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This volume is dedicated to Professor
Sreenivasa Rao Jammalamadaka.

Sreenivas has advanced statistical science

in deep and original ways, and the statistics
community has been enriched by his research
contributions, intellectual leadership, and
gracious mentorship of many

Jjunior colleagues.






Preface

Professor Sreenivasa Rao Jammalamadaka, formerly known as J.S. Rao and affec-
tionately known to most of us as JS, was born on December 7, 1944, at Munipalle,
India. Being under-aged for engineering studies was a blessing in disguise, and he
was among the first batch of students selected for the Bachelor of Statistics (B. Stat.)
degree at the Indian Statistical Institute (ISI), Kolkata. He received his Masters and
Ph.D. degrees also from the ISI, and has the distinction of being the first B.Stat.-
M.Stat.-Ph.D. of the ISI. He received his education from such legendary figures as
Professors P.C. Mahalanobis, J.B.S. Haldane, C.R. Rao, and D. Basu among others,
and worked with Professor C.R. Rao for his Ph.D. (1969) on path-breaking research
in the then newly emerging area of directional data analysis.

JS moved to the USA in 1969 and was a faculty member at the Indiana University
and then at the University of Wisconsin, Madison, before he moved to the Univer-
sity of California, Santa Barbara (UCSB) in 1976, where he has remained since
then. At UCSB he played a pivotal and leading role in creating the new Depart-
ment of Statistics and Applied Probability and was its first Chairman. He has been
a prodigious mentor to the graduate students in that department, having provided
guidance to as many of 35 Ph.D. students, at the last count. Throughout his career,
JS has been extraordinarily generous to his colleagues in India, inviting them to the
US and spending many of his sabbaticals helping Universities in India as well as in
other countries.

JS has published extensively in leading international journals. His research work
spans a wide spectrum which includes: goodness-of-fit tests, linear models, non-
parametric and semi-parametric inference, Bayesian analysis, reliability, spacings
statistics, and most notably, directional data analysis. He has written several books,
both for undergraduate students as well as for advanced researchers. He has collab-
orated with a large number of researchers from around the world in general, and
from India in particular. A Fellow of both the ASA and the IMS among other pro-
fessional organizations, he has served nobly the cause of statistics at many national
and international levels, including that of the President of the International Indian
Statistical Association.

The present volume consists of papers written by students, colleagues and collab-
orators of JS from various countries, and covers a variety of research topics which JS
enjoys and contributed immensely to. We are deeply thankful to these distinguished

vii



viii Preface

authors for their contributions. We thank the anonymous referees for their reviews
and suggestions. We also profoundly thank Dr. Kaushik Ghosh; his technical savvy,
editorial contributions, and deep dedication to this project was extremely impor-
tant in bringing this volume to fruition. Finally, it is a pleasure to acknowledge the
understanding, patience and cooperation of Springer-Verlag in bringing out this vol-
ume. It is with great pleasure and pride that we present to our readers this volume,
a Festschrift in celebration of the 65th birth anniversary of JS. We also take this
opportunity to wish JS many, many more years of active research which will enrich
the statistical sciences as well as foster further academic collaborations from far and
wide.

Ithaca, NY Martin T. Wells
Kolkata, India Ashis SenGupta
June 2010
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Chapter 1
Models for Axial Data

Barry C. Arnold and Ashis SenGupta

Abstract A variety of models have been proposed to accommodate data involv-
ing directional vectors in two-dimensions, with no identified start or end point. By
convention such directions are represented by points in the interval (0, ). Data
of this kind is called axial data. A survey of symmetric and asymmetric models
for axial data is provided here. In addition, for certain models, parametric inference
issues are addressed. In some cases, bivariate and multivariate extensions are readily
envisioned.

1.1 Introduction

To model directions of vectors which do not have an identifiable start point or end
point, an angular distribution with support (0, ) is required. It is customary that
such distributions are called axial distributions. Such axial data has often been ana-
lyzed by multiplying the observation values by two and then seeking a circular
distribution (often a circular normal density) that provides an acceptable fit. This
will be referred to as the “doubling” strategy, which is justifiable in many cases.
Some times alternative approaches can yield better results. An alternative approach
involves the recognition that in many cases axial variables can be viewed as circular
variables modulo 7. Arnold and SenGupta [1] proposed use of this approach, begin-
ning with a circular normal density for the original directions. Wrapping strategies
can be used to generate axial models as can approaches involving projections of
points determined by a given bivariate distribution in R2. In the case of circular data,
the most popular model is the von Mises or circular normal density. Its popularity
can perhaps be traced to the fact that the model is a two parameter exponential fam-
ily with minimal sufficient statistics that are simply sums of sines and cosines of the
observed values. Only by a doubling approach can a similar convenient exponential

B.C. Arnold (X)
Department of Statistics, University of California, Riverside, CA 92521, USA
e-mail: barry.arnold @ucr.edu

M.T. Wells and A. SenGupta (eds.), Advances in Directional and Linear Statistics, 1
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2 B.C. Arnold and A. SenGupta

family model be arrived at for axial data. This means that estimation and inference
for almost all axial models will be “computer intensive” to some degree.
It is time to begin the tour of “the world of axial modeling”.

1.2 Some Well-Known Circular Models

It will be useful to quickly review some of the more popular circular models (see
e.g., [6]). The first in our list (and undoubtedly the most frequently encountered) is
the circular normal distribution. A random variable ® will be said to have circular
normal distribution with parameters i € [0,27) and k¥ € (0, 00) if its density is of
the form

Jo(O; p. k) ocexple (6 — w)]I(0 < 6 < 2m). (1.1)

The parameter p corresponds to the mode of the density while « is a precision
parameter. If a random variable has density (1.1), we write &® ~ CN(u,k). An
alternative parameterization is possible, as follows

fo(8;a,B) xexplacosf + Bsin0]1(0 <6 < 2m). (1.2)

In this version the parameters o and B can take on any real value. Representation
(1.2), makes it evident that we are dealing with a two parameter exponential family
of densities and that, if we have a sample of size n from this density, the complete
minimal sufficient statistic will be

(Z:;l cos O, 27:1 sin @,-) . (1.3)

An alternative approach to the construction of models for circular data involves
“wrapping” a real variable or equivalently evaluating that variable modulo 27 to
yield a variable with support [0, 277). Thus we begin with a random variable X with
support (—oo, o0) and define the corresponding “wrapped” version of X by

® = X (mod 27). (1.4)

This is clearly a rich source of models since the distribution of X can be quite
arbitrary.

A popular choice for the distribution of the random variable to be “wrapped” is
a symmetric stable distribution. In particular, the normal distribution or the Cauchy
distribution is often selected. The wrapped normal distribution has a density express-
ible in the form of an infinite series, which complicates estimation and inference for
the model. On the other hand, a wrapped Cauchy distribution has a density of the
following relatively simple analytic form

fo@;a,B) o< (1 +acosf + Bsinf) 110 <6 < 2m) (1.5)
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where «, B are real valued parameters satisfying «? + 2 < 1. Note that the choice
a = 8 = 0 yields the uniform distribution on [0, 277).

Another distribution which is sometimes considered for univariate directional
data is the cardioid distribution with density of the form

fo(0;a,B) [l +acosf + BsinO]I(0 <6 < 2x) (1.6)

in which «, 8 are real valued parameters satisfying «?> + B2 < 1. A more general
model, introduced by [7] will be called a generalized Cardioid density. It is of the
form

fo@;a,B) <[l +acosf + Bsinf]I(0 <6 < 2x) (1.7)

in which «, 8, y are real valued parameters satisfying «? + 82 < 1. The third param-
eter y results in a flexible family of models which includes the wrapped Cauchy, the
uniform and the cardioid distributions as special cases correspondingto y = —1,0
and 1 respectively.

An alternative flexible family of circular distributions are those of the “angular”
form. For these , one begins with a arbitrary two-dimensional random vector (X, Y)
with support in the plane. We then consider the angle in the interval (0, 277) deter-
mined by the line joining the origin to the point (X,Y), ie.,, ® = tan"'(Y/X).
A popular choice for the distribution of (X, Y) in this construction is the classical
bivariate normal distribution.

The last entry in our list of general methods for creating flexible circular models,
is the technique of “restriction to the unit circle”. In this approach one begins with
an arbitrary joint density fo(x, y) whose support includes an open set that includes
the unit circle. Then one considers the values taken on by this joint density at all
points on the unit circle. Parameterizing the unit circle by 8 € [0, 27), one obtains
in this way a circular density of the form:

f(0) < fo(cosB,sinB)I(0 <6 < 2m). (1.8)

In fact, in this construction fy can be replaced by a quite arbitrary function of
two variables, not necessarily a joint density.

1.3 Introducing Asymmetry

Most of the circular models described in Sect. 1.2 are symmetric about their modes.
But of course, many real world data configurations do not exhibit such symmetry. To
accommodate this situation a variety of asymmetric versions of the densities have
been proposed. We will return to discuss asymmetry in more detail in the context
of axial distributions in Sect. 1.5. In the circular context, only a few brief comments
will be presented.
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First note that some of the constructions introduced in Sect. 1.2 clearly do not
always produce symmetric models. Thus, restriction to the unit circle and angular
constructions can easily yield asymmetric models. Likewise, wrapping an asym-
metric density usually leads to asymmetry (examples in the literature include the
wrapped exponential [5] and the wrapped skew-normal [9]).

Umbach and Jammalamadaka [10], motivated by [4] construction of skewed
distributions on the real line, describe a general method of constructing skewed
circular distributions. They begin with two symmetric circular densities f and g on
[—7, 7). Denote the distribution function of g by G and choose w to be an odd func-
tion with period 27 and satisfying |w(6)| < m. They then verify that the function
2 f(0)G(w(0)) is an asymmetric circular density.

Efforts to develop asymmetric versions of the circular normal distribution often
involve introduction of trigonometric functions of different frequencies. Thus, for
example, we might consider a model of the form

fo(0;a,B,y,6) xexplacosf + Bsinf + ycos(260) + §sin(30)]1(0 < 6 < 2x).
(1.9)

Analogous adjustments can be made to the generalized cardioid density to obtain
asymmetric versions.

1.4 Axial Models

As mentioned in the introduction, axial distributions are appropriate for modeling
the directions of vectors without an identifiable beginning or end point. They typ-
ically will take on values between 0 and . Two general strategies are considered
for generating axial models from available circular models: wrapping and doubling.
The doubling approach involves multiplying the axial variable by 2 and assuming
that the doubled variable has one of the available circular distributions described in
Sect. 1.2. Thus if our axial random variable is denoted by ® and if g(0) is a given
circular density, then the density for ® will be of the form

fo(0) =2g(20)1(0 <0 < ). (1.10)

Generally speaking such models will be suspect, because there seems to be little
reason to expect that two times an axial variable will be sensibly modeled by a well
known circular distribution. Nevertheless this technique does provide a very wide
ranging flexible array of possible models which might be useful for modeling and/or
prediction. There is an exceptional case in which doubling does seem to make sense,
to be described later.

The second approach for generating axial models involves wrapping, but in this
case wrapping around a half circle instead of a full circle as was done to generate
circular models. Thus one begins with an arbitrary real valued random variable X
and defines an axial variable ® by setting



1 Models for Axial Data 5

® = X (mod ). (1.11)

Generally speaking it is desirable that axial density f(0) should have the property
that limg_,, f(6) = f(0), since the angles 0 and 7 are indistinguishable. In parallel
fashion it is reasonable to expect that a circular density will satisfy limg_,,, f(6) =
£(0). Such restrictions will limit the class of acceptable densities for the random
variable X that is to be wrapped.

Perhaps the most natural genesis for an axial variable involves envisioning a
circular random variable that is imperfectly visible in the sense that any pair of
values of the form 6 and 8 + 7 are indistinguishable. Thus if the original circular
variable is denote by @ the density of the observed axial variable will be of the form

Jo(0) =[fe(0) + fo (0 + )] 1(0 <0 < 7). (1.12)

An alternative viewpoint, is that this density results from the fact that ® =
@ (mod ).

A particularly popular version of this construction begins with circular normal
distribution for @ and results in what is called an axial normal distribution for ® of
the form

fo(0) «x cosh(asinf 4+ BcosO) I(0 <6 < ). (1.13)

See [1] for more detailed discussion of such axial normal variables. These axial
normal variables, derived in a natural fashion from circular normal variables, are
somewhat more difficult to deal with because the corresponding collection of
densities does not comprise an exponential family.

In passing, it may be noted that beginning with a circular normal density and
using the doubling approach does lead to an exponential family of axial densities of
the form

fo(0;a, B) x explacos20 + Bsin20] I(0 < 6 < m). (1.14)

The attraction of such models, with their conveniently available minimal sufficient
statistic, is undeniable. Despite the lack of logical justification for use of the dou-
bling approach in this context, it must be admitted that they comprise a flexible two
parameter family of axial distributions which merits consideration for use in fitting
data.

Instead of using the wrapping or the doubling approach, angular axial models
may be considered. To this end we, as in the circular case, begin by considering a
bivariate random vector (X, Y) with support in the plane. Then let ® denote the
angle in the interval (0, ) determined by the extended line joining the origin to
the point (X,Y). Thus @ = tan~!'(Y/X) + (r/2) where tan™! is chosen to range
over the interval (—m/2,7/2) as is customary. Although the model is motivated
by consideration of angles determined by a random point in the plane, it can be
described in a slightly simpler fashion. The random vector (X, Y) only enters into
the model in terms of its ratio ¥/ X and since the distribution of (X, Y) is arbitrary,
so is the distribution of the real random variable W = Y/ X. So the general angular
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axial model can be represented in the form
O =tan ' W + (/2) (1.15)

where W is an arbitrary real random variable.

A popular choice for the distribution of (X, Y) in this construction is the classical
bivariate normal distribution with mean vector i and variance covariance matrix X'
With this choice of distribution for (X, Y) the random variable W = ¥/ X will have
a Cauchy distribution and as u and X' range over all possible values, the location
and scale parameters of the Cauchy random variable W will range over all possible
values, i.e., —00 < i < oo and 0 < 0 < oo.

With such a choice for the distribution of (X,Y), or equivalently of W, the
resulting axial distribution is known as the angular central Gaussian distribution [8].
There are several possible parameterizations but the one providing the most simply
interpretable parameters is of the form

fo() o (1+asin (z (9+%—ﬂ)))_1 10<6<m).  (1.16)

The parameter o controls the precision (or peakedness) of the density while S
controls the location of the mode.

One obvious advantage of this model is that a simple transformation applied to
the density yields a Cauchy density so that inference techniques appropriate for
Cauchy distributed data may be directly used in analysis. But there is another,
perhaps unexpected special property of the angular central Gaussian (ACG) dis-
tribution. These features of the model become more apparent when alternative
parameterizations are considered.

A parameterization of the ACG density that highlights its linkage with a
Cauchy(u, o) density is as follows:

20 I(0<0 <)

. 2 —g2
7(l+0%+ pu?)[l - 1+022—M+u2 sin(26 — ) — ;-FZT-F(:LZ cos(26 — m)]
(1.17)

Jo(0) =

where —oo < 4 < ocand 0 < 0 < 0.
Instead one might consider a version of the ACG density parameterized as

follows
V1=82—y2 I(0<0 <m)

Jo(0) = (1 + ysin(20) + § cos(26))

(1.18)

where the parameters y and § are constrained by
0<y>+6%<1.

Of course the choice y = § = 0 leads to a uniform distribution on [0, 7). If ® has
the density (1.18), it is clear that the doubled random variable A has a density of
the form
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Fay(d) o< (14 ysind +8cos )™ 1(0 < A < 27), (1.19)

which is recognizable as a wrapped Cauchy distribution (c.f., (1.5)). It would thus
be possible to double data arising from an ACG distribution and analyze the doubled
data using a wrapped Cauchy model.

Remark. The double linkage between the ACG distribution and the Cauchy distri-
bution raises an interesting characterization problem. It is apparent from the above
discussion that if W has a Cauchy(0, 1) distribution, then for some a, b the random
variables U = a +bW (mod 27r) and V = 2(tan" ! W + (n/2)) = 2tan ' W +
are identically distributed. Is this only true in the case in which W has a Cauchy
distribution?

Parallel to the development of the generalized cardioid circular model, it is natu-
ral to extend the ACG distribution by introducing an additional parameter, say p, to
consider generalized ACG densities of the form

fo(0) < [1 + ysin(20) + §cos(26)]° 1(0 <0 < 7). (1.20)

Yet another manner in which an axial density can be constructed is by “restriction
to the unit half-circle”. As in the analogous method of generating a circular density,
one begins with an arbitrary joint density fo(x, y) whose support includes an open
set that includes the unit half circle. Then one considers the values taken on by this
joint density at all points on the unit half circle. Parameterizing the unit half circle
by 8 € [0, ), one obtains in this way an axial density of the form:

f(0) x fo(cosB,sinB)I(0 <6 < ). (1.21)

1.5 Asymmetric Axial Models

For axial distributions, as was the case for circular models, the majority of the
popular densities are symmetric about their modal value. Asymmetric variations
are however available. Wrapping an asymmetric real random variable X around
the unit half circle will typically yield an asymmetric axial distribution. Such is
also the case if we make use of the angular projection approach, i.e., consider
©® = tan~!' W + (/2) where W is an arbitrary real random variable. In this con-
struction, the choice of a Cauchy distribution for W will lead to a symmetric density
for @, but other choices for the distribution of W lead to asymmetric axial densi-
ties. Even a standard normal choice for W leads to asymmetry! An intriguing open
problem is to characterize the class of distributions for W that will lead to symmet-
ric axial distributions. It is not difficult to verify that this class includes t-densities
with possibly fractional degrees of freedom, but presumably there are many more
possibilities.

Finite mixtures of symmetric axial densities will typically exhibit asymme-
try, as will finite logarithmic mixtures. More generally, if f1(0), f2(8),..., fm(6)
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are m symmetric axial densities, then subject to non-negativity and integrability
constraints one may consider the following usually asymmetric axial models

f(6) [Z a; f; (9)} JI(0<6 <), (1.22)

i=1
and
m
1) « [H{ﬁ- (9)}“1} /100 <6 <). (1.23)
i=1
It is possible to develop asymmetric versions of the axial normal distribution

by the introduction of trigonometric functions of different frequencies. Thus, for
example, one might consider a density of the form

fo(8;a,B,y,8) « cosh[asinf + B cosO + ysin(20) + § cos(30)]1(0 < 0 < )
(1.24)

Analogous adjustments can be made to the ACG and extended ACG model to obtain
asymmetric versions such as

fo(0) < [1 + ysin(20) + 8 cos(26)) + A sin(40) + t cos(66)]° 1(0 < 0 < 7).
(1.25)

1.6 Bivariate Axial Distributions

Bivariate (and multivariate) versions of the axial distribution (1.15) are readily
constructed by choosing (W;, W5) to have a bivariate (or W to have a multi-
variate) Cauchy distribution with Cauchy(0, 1) marginals and then applying a
transformation of the form (1.15) to each coordinate, i.e., define

O; = tan ' (uy + o1 Wy) + (r/2)

for eachi.
For some applications it may be desirable to have a bivariate (or multivariate)
axial distribution with conditional densities belonging to specific axial families.
For example, if we wish to deal with bivariate distributions with conditionals in
the generalized ACG family (1.20), we may consider densities of the form

fo,,0,(01,62) x [(1, sin 2601, cos 2601) A(1, sin 26;, cos 292)’]p
1(0<6; <7 0<6,,m). (1.26)
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Itis evident that (1.1) does indeed have all conditional densities of ®; given ®, and
of ®; given O in the family (1.20). The choice p = —1, yields densities with ACG
conditionals.

Similarly, it is possible to describe a bivariate density with all conditionals in the
axial normal family (1.13). It will be of the form;

f(61,65) o cosh[(sin 1, cos 01)B(sin 85, c0s 62))1 1(0 < 0; < 7,0 < 6, < ),
(1.27)
where B is a 2 x 2 matrix of parameters.
In the case of axial model (1.14), the fact that this is two parameter exponential
family allows us (using [3]) to identify the class of all bivariate axial densities with
conditional densities of the form (1.14). Such densities will be of the form

f(61,62) = exp][(1,sin 201, cos 261)A(1, sin 265, cos 26,)']
1(0< 6y <m,0<6,,m), (1.28)

where A is a 3 x 3 real matrix in which a;; is a normalizing constant that is a
function of the other a;;’s so that the density will integrate to 1. Multivariate versions
of this construction can be analogously defined.

Asymmetric multivariate axial models can be constructed using basically the
same devices that were used to construct asymmetric univariate axial models. More
details on these constructions may be found in [2].
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Chapter 2

Asymptotic Behavior of the Universally
Consistent Conditional U-Statistics

for Nonstationary and Absolutely Regular
Processes

Echarif Elharfaoui, Michel Harel, and Madan L. Puri

Abstract A general class of conditional U-statistics was introduced by W. Stute
as a generalization of the Nadaraya—Watson estimates of a regression function. It
was shown that such statistics are universally consistent. Also, universal consis-
tencies of the window and kj,-nearest neighbor estimators (as two special cases
of the conditional U-statistics) were proved. Later, (Harel and Puri, Ann Inst Stat
Math 56(4):819-832, 2004) extended his results from the i.i.d. case to the absolute
regular case. In this paper, we extend these results from the stationary case to the
nonstationary case.

2.1 Introduction

Let {Z; = (X;,Y;); i € N*} be a sequence of random vectors with continuous
distribution functions H;(z), i € N*, z € R? x R®, defined on some probability
space (£2, A, P).

Assume that H; admits a strictly positive density and H; has the two marginals
F; and G;.

Let /1 be a function of k-variates (the U kernel) such that forsomer > 2,h € L},
which means that E{supg |h(Yp)|"} < +o0 (where sup extends over all permuta-
tions B = (B1,...,PBr) of length k, that is, over all pairwise distinct f1, ..., Bx

taken from N*) which implies that for all integers i1, i2,...,if (i1 < iz < ... <
ix) h(Y;,,..., Y ) € Ly, the space of all random variables Z for which |Z|" is
integrable. In order to measure the impact of a few X’s, say (Xy,..., Xg), on a
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function A(Y1, ..., Yy) of the pertaining Y’s, set
m(x) = m(xy,...,xx) := E[h(Y1,...,Y)| X1 = x1,..., X = x¢] 2.1

where m is defined on R%¥.
For estimation of m(x), [7] proposed a statistic of the form

Yph (¥, ... . Yg) TT5—y KI(x; — Xp,)/ hnl
Y g [T52 Kl(xj — Xp,)/ ha]

un(X) = up(x1,...,X5) = 2.2)

where uy, is defined on R%* K is the so-called smoothing kernel satisfying [ K (u)
du = 1 and {h,, n > 1} is a sequence of bandwidth tending to zero at appropriate
rates. Here summation extends over all permutations 8 = (81, ..., Br) of length k,
that is, over all pairwise distinct 81, ..., B taken from 1, ..., n. Stute [7] proved the
asymptotic normality and weak and strong consistency of u, (x) when the random
variables {(X;, Y;), i > 1} are independent and identically distributed. Harel and
Puri [3] extended the results of [7] from independent case to the case when the
underlying random variables are absolutely regular. Stute [9] also derived the L,
convergence of the conditional U -statistics under the i.i.d. set up.

If a number of the X;’s in the random sample are exactly equal to x which can
happen if X is a discrete random variable, PY (-|X = x) can be estimated by the
empirical distribution of the Y;’s corresponding to X;’s equal to x. If few or none
of the X;’s are exactly equal to x, it is necessary to use Y;’s corresponding to X;’s
near x. This leads to estimators ﬁ,{ (-|X = x) of the form

n
PYCIX =x) =) Wai(0)lly, e

i=1

where W, (x) = Wyi(x, X1,...,X,) (1 < i < n) weights those values of i for
which X; is close to x more heavily than these values of i for which X; is far from x
and 114 denotes the indicator function of A.

Let g be a Borel function on R* such that g(Y) € L,. Corresponding to W, is
the estimator /,, (x) of [(x) = E(g(Y)|X = x) defined by

In(x) = Z Wai(x)g(Yi).

i=1

More generally if we now consider the estimates of m(x) defined in (2.4), this leads
to weighting those values of 8 for which Xg = (Xg,,..., Xp, ) is close to x more
heavily than the values of 8 for which Xg is far from x.

This is why, as in [8], we study a fairly general class of conditional U -statistics
of the form
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ma(x) =Y Wpu(x)h(Yp) (2.3)
B

designed to estimate m(x), where Wg ,(x) is defined from a function W, (x,y) by
Wgn(x) = Wu(x,Xg), Yg = (Yp,,...,Yg,), and the summation in (2.3) takes
place over all permutations 8 = (B4,...,Px) of length k such that 1 < ; <
n,i=1,...,k.

Remark 2.1. The estimator defined in (2.2) is a special case of the estimator defined
in (2.3), see (2.23).

In order to make m, (x) a local average, Wpg , (X) has to give larger weights to those
h(Ypg) is close to x. For this general class of conditional U -statistics (defined in
(2.3)) and for i.i.d. random variables, [8] derived the universal consistency. Harel
and Puri [4] extended his results from the i.i.d. case to the absolute regular case.
In this paper, we extend it to the nonstationary case and absolutely regular r.v.’s
which allow broader applications that include, among others, hidden Markov models
(HMM) described in detail in [4].
We shall call Wg ,, universally consistent if and only if

my(X) > m(X) in L,

under no conditions on % (up to integrability) or the distribution of {(X;, ¥;), i > 1}.
Here X=(X?,..., X,?) is a vector of X ’s with the same distribution as (X1, ..., X%)
and independent of {(X;, Y;), i > 1}.

2.2 Preliminaries

Let (Z;);>1 be a stochastic process indexed by the positive integers, taking value
in a finite dimensional Euclidean space H. Identifying H with a product of a finite
number copies or the real line, we write H; for the distribution function of Z;. We
will assume that the process has some form of asymptotic stationarity, implying that
the sequence H; converges in a sense to be made precise to a limiting distribution
function H. _

Fori < j, let .A{ denote the o-algebra of events generated by Z;, ..., Z;. We
shall say that the nonstationary stochastic process is absolutely regular if

sup max E { sup |P(A|.A{)—P(A)| =Bk)* L0 as n — o0

1<j<n—k
neN* 1=j=n Ae.A‘j?°+k

where N* = {1,2,...}.
All along the paper, we assume that (*) holds with a geometrical rate;
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Z mﬂlsﬁ(m) < oo forsomed > 0. (2.4)

m>1

We consider a parameter £ in H whose components can be naturally estimated by
U-statistics. To be more formal and precise, we assume that £ is defined as follows.
Let k be an integer, to be the degree of the U-statistics. Let ¢ be a function from HF
into HI, invariant by permutation of its arguments. We are interested in parameters
of the form

k
= dH®* = SO dH(z). 2.5
e=[ o IRE w [Jart @5

and the function ¢ is called the kernel of the parameter &.

Example 2.1. Take H to be R. The mean vector corresponds to taking k = 1 and ¢
is the identity.

Example 2.2. Take H to be R2. Consider £ to be the two-dimensional vector whose
components are the marginal variances. We take k = 2 and ¢ is going to be
a function defined on (Rz)z. It has two arguments, each being in R2, and it is
defined by

2 2

u? +u' L VY ,

, -w].
2 2

¢ ((Lt, V)’ (ulv V/)) = ( — uu

Such a parameter can be estimated naturally by a U-statistics, essentially replac-
ing H®* in (5) by an empirical counterpart. By using the invariance of ¢, the
estimator of £ is then of the form

-1
£, = (Z) > $(Zp,..... Zg)- 2.6)
p

To specify our assumption on the process, it is convenient to introduce copies
of H. Hence we write H;, i > 1, an infinite sequence of copies of H. The basic idea
is to think of the process at time i as taking value in H; and we think of each Hj; as
the ith component of H*. We then agree on the following definition.

Definition 2.1. A canonical p-subspace of H* is any subspace of the form H;, &
... ®H;, with 1 <iy <--- <i,. We write S, for a generic canonical p-subspace.

Remark 2.2. For (iy,....ip) # (j1.....jp) if wenote S, = H;, &... ®H;, and
S, =H;, &... H;,, wehave S, # S/, with S, C H* and S, C H*.

The origin of this terminology is that when H is the real line, then a canonical
p-subspace is a subspace spanned by exactly p distinct vectors of the canonical
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basis of H. We write } 5 for a sum over all canonical p-subspaces included
in H".

To such a canonical subspace S, = H;, & ... H; , We can associate the distri-
bution function Hs, of Ziy,.... Z; p) as well as the distribution function with the
same marginals

H®? = ®i<j<pH;, = Qu,cs, Hi. 2.7)

i

Clearly the marginal of H ®S? are independent, while that of Hs , are not.

Consider two nested canonical subspace S, and Sg_, where Sx_, C H" ©S,,.
For a function ¢ symmetric in its argument and defined on S, &Sk _ ,, we can define
its projection onto the functions defined on S, by

L€S) = (2 Sk_p) = /S B, y) AHDS1 (). 2.8)

Identifying S, ® Sk—, with H* and H? with S p» that allows to project functions
defined on H¥ onto functions on HPZ. However, with this identification, the projec-
tion depends on the particular choice of Sg_, in H". To remove the dependence in
Sk—p, we sum over all choices of Sy, in H" © S by

-1

n—

s, (2) = (k _ p) Yo @Sy (2.9)
p Sk—pCH'OS)

Let k be an integer and for each n > k, consider a kernel ¢, = ¢ of degree k

depending on n.
A U-statistics of degree k is defined by

-1
Uy = (Z) > $a(Zpyo . Zp)s (2.10)
5

we can then define an analogue of Hoeffding decomposition when the random vari-
ables come from a nonstationary process. For this purpose, consider, firstly, an
expectation of U, if the process had no dependence, namely,

-1
Upo = (Z) S | ¢ dH®x. @2.11)
Sy CH”? Sk
Then forany p = 1,..., k, we define
-1
n
Un,p = ( ) > [ $s, d ®s,cs, 6z, — Hy) (2.12)
PJ) s, cun /e

where 4y ; is the Dirac function.
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Finally, for p > k, we set
Un,p =0. (2.13)

The analogue of Hoeffding decomposition is the equality

Un= Y <k>U,,,,,. (2.14)

0<p<k P

Note that this decomposition makes an explicit use of convention (2.13), and this is
why this convention was introduced.

We now need to specify exactly what we mean by asymptotic stationary of a pro-
cess. For this, recall the following notion of distance between probability measures.

Definition 2.2. The distance in total variation between two probability measures
P and Q defined on the same o -algebra A is

|P — Qla = sup |P(4) — Q(A4)].
AcA

If S, is a canonical subspace of H*, we write o5, the o-algebra generated by the
Z;’s with H; C S,. We write P the probability measure pertaining to the process
(Z;)i>1, which is a probability measure on H*.

Definition 2.3. The process (Z;);>1 with probability measure P on H> is geo-
metrically asymptotically stationary if there exists a strictly stationary process with
distribution Q on H®°, and a positive t less than 1, such that fori > 1,

|P = Qloy, <7 (2.15)

We suppose that there exists a strictly stationary process (Z);>1 with probabil-
ity measure Q on H, which is absolutely regular with the same rate as the process
(Zi)i=1. H is the distribution function of Z, H admits a strictly positive density
and H has the two marginals F and G.

We define the function ¢* on H; by

zeH; — ¢*(z HF 0 H,)) = / ¢z, y) dH®*D, (2.16)
HK OH;

Next, we denote

n
Ur =n" Z/H ¢*d(zx — H).
1

i=1

2.3 Assumptions and Main Results

In this section, we identify H = H’' x H” with R? x R. For a generic canonical
p-subspace S, of H*®, we write S; x and S, x its projections respectively in H'*®
and H"*°.



2 Asymptotic Behavior of the Universally Consistent Conditional U-Statistics 17

We consider the nonstationary sequence of random vectors {Z; = (X;,Y;); i €
N*} with values in R4 x RS and continuous distribution functions H; and H; has
the two marginals F; and G;.

We assume that the sequence {Z;};>1 is absolute regular with rates (2.4) and
(2.15) is satisfied with its associated stationary sequence {Z = (X Y ); i € N*}
of stationary random vectors.

For the ease of convenience, we shall write Wg for Wpg ;.

Consider the following set of assumptions:

(i) There exists functions Vj(x,y) on R24¥ such that for each [ € Lcr, 2™ =
(2152 20) € R and y™ = (y1,..., yy) € R*"

2.p Vn(x.2p)l(yp)
> p Va(x,2p)

Z Wy (X’ Zﬁ)l(yﬂ) =
B

where zg = (z8,,...,28,) and yg = (¥g,, .., Vg )-
(ii) There exists a function V(x) on R4¥ such that for each scalar function q on R4k
verifying

/WﬁM@WﬂﬁW@<m

we have

-1
lim (Z) Z [ q(2)Vy(x,2)dF®S1% (z) = q(x)f(x)/ V(z)dz

n—o0
S1.xCH"

where f (x) = ]_[];-:1 f(x;)and f is the density function of F.
(iii) Define the kernel of degree k by

$r(23) =B Va(x.2) | [ Valx P ),

Suppose that
sup [ |pul*T? dPoy, < 00 2.17)
Sk CH®® JSg
sup [ |pa*T dQy, < o0 (2.18)
Sk CH®® JSg
where § > 0.

Remark 2.3. Our conditions (i) and (ii) are completely different from conditions
(ii) to (v) in [8]. Our conditions are more general and more easy to verify. More, the
condition (i) in [8] is not necessary.

The following theorems generalize Theorems 2.1, 2.2, 2.3 and 2.4 in [4] from the
stationary dependent case to the nonstationary dependent case.
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Theorem 2.1. Assume that h € L. Then under (i)—(iii), (2.4) and (2.15),
my(X) > m(X) inL,, asn — o0,
where r = 2 + 26, that is
E [|my(x) — m(x)|" u(dx)] > 0asn — oo (2.19)

where 1 denotes the distribution of (X1, X2, ..., Xg).

Corollary 2.1. Under the conditions of Theorem 2.1 and (2.34) in Sect. 2.4, my(X)
— m(x) with probability one for p-almost all x.

Remark 2.4. In [4], we supposed that & is bounded, this condition is not necessary
now.

Theorems 2.2 and 2.3 deal with two special cases: window weights and NN-weights.
Consistency of window estimates for the regression function has been obtained
by [2] and [5]. NN-weights for the regression function have been studied in [6],
Theorem 2.

In what follows, | - | denotes the maximum norm on R¢. We also write

Xs — x|| := max |Xg — x;|.
IXp — x| = max |Xp, —xil

To define window weights, put (see [8])

Wp(x) = (1)1[||Xﬁ—x||shn]/ 2_p Miixg—xl<hs)  if well defined (2.20)

otherwise.

Here h, > 0 is a given window size to be chosen by the statistician. Then we have
the following results:

Theorem 2.2. Assume h,, — 0 and nh,“l’ — oo as n — oo. Then, under the
conditions (2.4) and (2.15), we have

my(X) > m(X) in L,,

where Wg(X) in (2.3) is given by (2.20).

For the NN-weights, recall that X ; is among the k,NN of x € R? if d;(x) :=
| X; — x| is among the k,-smallest ordered values d.,(x) < ... < du:n(x) of the
d’s. Ties may be broken by randomization.

For a given 1 <k, <n, set

k;d if Xg. is amongthe k, — NN of x; for1 <i <k

W, =
p(x) 0 otherwise.

2.21)
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Theorem 2.3. Assume that k, — oo and k,/n — 0 as n — co. Then, under the
conditions (2.4) and (2.15), we have

my(X) > m(X) in L,
where Wg(X) in (2.3) is given by (2.21).
We now consider as estimator of m(x), the statistics of the form
My (X) = utn(X) (2.22)

where u, (x) is defined in (2.2). Then, in view of (2.3) we have

151 Kl(xj — Xp,)/ ha]
> TT5—1 Kl(x; — Xp,)/ ]

Wp . (X) = (2.23)

where K(x) is a so-called smoothing kernel satisfying [ K(u)du = 1 and
limy—oo [u|K(u) = 0 and {h,, n > 1} is a sequence of bandwidths tending to
zero. This special case was studied by [7] for i.i.d. random variables, and from
Theorem 2.1, we can generalize his result for nonstationary dependent random vari-
ables. The following theorem establishes that the universal consistency still holds
for conditional U-statistics involving kernel K and a sequence of bandwidth /,,.

Theorem 2.4. Assume that h,, — 0 and nhg — oo asn — o0o. Then, under the
conditions (2.4) and (2.15), we have

my(X) > m(X) in L,,

where m(x) is given (2.1).

2.4 Proof of Theorems and Corollary 2.1

First, we show that m,, is the ratio of two U -statistics. Let x = (x1, ..., x¢) be fixed
throughout. Let

-1
Un(h,x) = Up(x) = U, = (Z) > h(Yp)Vax, X,g)// Vo (x, u)dF®* (u).
5

Hence m, (x) = Uy (h, X)/U,, (1,x) and U, (h, x), for each n > k, is a nonstationary
U -statistic as defined in (2.10) with a hind depending on 7.
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Consider the sequence of functionals

—1
uhx)=0,= (") Y [ puar®%
k S, chn Y Sk
k

where ¢, is defined in (iii).
Note that 68, = E(U,).
The decomposition defined in (2.14) can be written as

“ (k
U”:9"+Z( )Un,p
=1 \7

where U, p is defined as in (2.12).
To prove Theorem 2.1, the following lemmas are needed.

Lemma 2.1. Under the conditions of Theorem 2.1
E(Un,p)* = O(n™?).
Proof. We shall consider the case p = 2. The proofs in the cases ¢ = 3,...,k

are analogous and so they are omitted.
We first note that

-1
n
Un,2 = (2> Z / ¢52d®]}-]1ics2 (82,- - Hl')
S, CH” S2
so we have
-1
n
E(Uyp)* = (2) Z Z J((i1,12), (I1,12))
1<iy<iz<mn 1<ly<lr<n

where
J((i1,i2), (I, 1)) = E (/S ¢S2d®15,-52H,-j (SZl.j —H;)
2
x/ bs;de| <oy, 8z, — Hlm))
S5
S> = H;, ® Hj, and S; = H;, & Hj,.

So from condition (2.4) and condition (iii), we have from Lemma 2.1 in [10] the
inequalities:
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DOIf1 <iy <ip <l <l then

J((r.i2). (. 1)) < M{B(la — 1)}T98: Iy — Iy > i — iy

and
J((i1.i2). (11, 12)) < M{B(ia — i)} T8 iy —iy > o — Iy

where M is a finite positive constant.
Thus, using (2.24) and (2.25), we obtain

> J((i1.i2). (. 1)) | = O(n?).

1<iy<ipz<li<lz<n

Similarly
i) If1 <iy <ly <ip <ly <n,then

> J((i1,i2), (I1,12))| = O?).
1<iy<ly<iz<lr<n
G If1 <iy <y <1, <ir <n,then
Yo JlGhi2), (k)| = O(?).
1<ii<li<lx<iz<n
From (2.26), (2.27) and (2.28), we obtain
E(Un2)? = 0(n™?).
Thus the result for the case p = 2 is proved.

Lemma 2.2. Under the condition of Theorem 2.1, for p-almost all x

On(h,x)
On(1,x)

— m(x), n — oo.
Proof. By definition, we have

n

-1
O (h,x) = 0 = (k) > PndH®SK
Sy CH" Sk
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(2.24)

(2.25)

(2.26)

(2.27)

(2.28)
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Put .
6! (h.x) = (Z) / h(y)Vy (x, 2)dF®5% (y, z).
S CH”
From condition (2.15), we deduce
|6, (h,x) — 6, (h,x)| > 0 asn — oco. (2.29)

From condition (ii) in Sect. 2.3, we have

Jim (Z) Z /S m(z) Vi (x, 2)dF®S1% (z) = m(x) f (x) / V(z)dz

K CH”

(2.30)

and so

Tim_ (Z) ZCJHI’” [S Vo (x, 2)dF®S15 (z) = f(x) / Vz)dz. (2.31)

By definition, we have

en<h,x)=<2) > h(y)vn<x,z)dF®Sk<y,z)

SCH
) (”)_ Z/s (/S” E(h(y)|X = 2)Va(x, Z)dG®S2,k(y)>
xdF®ka2H;
: <Z)_l z [ mEVa o). (2.32)
§1 4 CHI"

From (2.29)-(2.32), we deduce easily that

0n (R, x)
0,(1,%)

— m(x), n — oo.

To prove Theorem 2.1, from Lemmas 2.1 and 2.2, we now have to show that for
p-almost all x,
Uy.,1(h,x) — 0 in probability.
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Since
Unp(h,x) =n"" Z ¢s,d ®m;cs, 6z, — H;)
S| CH” S1
n
=n"! Z/ ¢H,‘d(82i — H;)
i=1"Hi
we have

n 2
W) =2 (87, — H;
EUn1)? = n E(Z[Hi"bﬂ' 5z H))

i=1

n 2
= -2 d 8 . i
n ;E([Hi@}ﬂ, 0z H))
+ 2n~2 Z E (/ ¢Hid(82i - Hl))
H;

1<i<j<n

x( ¢, d(8z; —Hj))} )
H

From Lemma 2.1 of [10] and condition (iii), we have

E(Un,1)* <2n7nM(2,h) + 4’1_2M‘7}F’S(V7h) Xn:(P + 1),3%(17)
p=1
—om™)
where M (t,h) = E{supg |¢(Xp. Yp)|'}, which implies
E(U,1)*=0m™). (2.33)

From Lemmas 2.1 and 2.2 and from (2.33), we have

Uy, (h,x) — m(x)f(x)[ V(z)dz

and
Un(1,x) > f(x) / V(z)dz in probability

as n — oo for w-almost all x.
It remains to prove the uniform integrability.
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It is an easy convergence of the Jensen’s inequality

r

sup E 1| > Va(X.Xp)h(Yp)l/ Y Va(X. Xp)
B B

neN*

< sup £ D Ve Xp)h(Yp)I"/ D Va(X. Xp)
neN* 8 8

< E {sup|h(Yp)|"p < +oc.
B

and Theorem 2.1 is proved.
The proof of Corollary 2.1 is a consequence of Lemma 2.1 and Lemma 2.3 below.
For a d-dimensional vector V', consider the norm || V|| = max;<;<g | V)|, This
norm is equivalent to the Euclidian norm and easy to work with here. We will use
this norm in Lemma 2.3 below see also [1].

Lemma 2.3. Let (V,)n>1 be a sequence of d-dimensional centered absolutely
regular and non necessarily stationary random vectors with rate satisfying

Y () B < oo (2:34)
i>1
suIIDE(IIViII’) < oo. (2.35)

Then .
n! Z Vi — 0 with probability 1, as n — oco.

i=1
)

> e) . (2.36)

) . (2.37)

Proof. For € > 0,

P(wl=e) = 7 (ms,
< Y P(

1<j=<d

n

1 .

i=1

1 (;

i=1

Forall 1 < j < d, one has from Markov’s inequality that

1
e'n’

n

EE:IGU)

i=1

o (;

i=1
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By Lemma 5.2 of [3], one has that

n r
E ( Pk ) <cn'l? (2.38)

i=1
From the above two inequalities, one deduces that

n

,
- C
ElD_ v ) < =nl2 (2.39)

=
i=1

Since r/2 > 1, the last inequality implies that forall 1 < j <d,

n
ZP(%ZVI-U) ze><oo

n>1 i=1

which, in turn, implies that

n
ZP %ZVI >e | < o0.

n>1 i=1

Lemma 4.3 then follows by Borel-Cantelli theorem.

The proofs Theorems 2.2 to 2.4 are also consequences of Theorem 2.1 by using
technics similar as in the proofs of Theorem 2.2 to Theorem 2.4 in [4]: that is to
verify that conditions (i)—(iii) are satisfied.
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Chapter 3

Regression Models with STARMA Errors:
An Application to the Study of Temperature
Variations in the Antarctic Peninsula

Xiaoqi Gao and T. Subba Rao

Abstract Motivated by spatio-temporal problems that occur in many areas such as
environment, geography etc., we propose multivariate regression models with space-
time ARMA errors. The methods of model identification and estimation are studied.
The asymptotic properties of the estimators have been derived and simulations are
provided. The methodology is applied to the analysis of monthly mean surface tem-
peratures at five locations in the Antarctic Peninsula. This area of Antarctic is of
great concern to climatologists, as it is believed that there is higher rate of warming
compared to the rest. During the period from January 1978 to December 1998, the
temperatures at all the five locations in the Antarctic Peninsula have increased. Sub-
stantial warming were detected at Faraday/Vernadsky and Rothera stations with the
warming rate of 1.07 °C/decade and 1.08 °C/decade respectively and both trends are
significant at level 1%.

3.1 Introduction

The Intergovernmental Panel on Climate Change (IPCC) confirmed that the global
average surface temperature has increased by 0.61+0.2 °C since the late 19th century
(see [4]). Though the earth is unequivocally warming, the geographical patchiness
of the recent climate change is evident since it has been reported that the Antarc-
tic Peninsula (the northern most part of the mainland of Antarctica) is one of the
rapid warming areas in recent 50 years (see [4]). Since the temperature records
at industrial areas may be masked by both the increased concentrations of short-
lived sulphate aerosols and the heating of urban meteorological stations, it has
been suggested by [13] that the temperature trends of Antarctic stations are a par-
ticularly important indicator of climate change. Furthermore, the warming of the

X. Gao (X))

Mathematics Department, University of Manchester, Alan Turing Building, Oxford Road,
Manchester, M13 9PL, UK

e-mail: Xiaoqi.Gao@postgrad.manchester.ac.uk

M.T. Wells and A. SenGupta (eds.), Advances in Directional and Linear Statistics, 27
DOI 10.1007/978-3-7908-2628-9_3, (© Springer-Verlag Berlin Heidelberg 2011


Xiaoqi.Gao@postgrad.manchester.ac.uk

28 X. Gao and T.S. Rao

coastal areas in the Antarctic will lead to a collapse of ice shelves which results
in an increase of sea level. Therefore, the analysis of temperatures at the Antarctic
Peninsula is critical for understanding the dynamic behavior of climate change.

There are a number of recent studies concerned with the temperature trends of
the Antarctic Peninsula (see, e.g., [12, 13]). These studies agree that the Antarctic
Peninsula is warming and the statistically significant warming happened at Fara-
day/Vernadsky and Orcadas stations with the rates of 0.5 °C/decade and 0.2°C/
decade respectively. These trends were obtained by fitting a simple least square
regression model with independent errors. Since the temperature series is obvi-
ously temporally autocorrelated, the significance of the trend using the adjusted
t-test [9, 11] is not appropriate.

It is widely believed that besides temporal autocorrelation in the temperatures
at each location, there is a strong correlation between spatial locations (see [5]). In
time series modeling, these facts need to be taken into account. In view of this, the
temperatures at different locations in the Antarctic Peninsula is obviously a spatio-
temporal problem. Based on the belief that we will have a better insight into the
dynamics by including spatial information in modeling rather than analyzing each
series separately, we propose a multivariate regression model with correlated errors
having spatio-temporal structure which takes into account both spatial dependen-
cies and temporal autocorrelations. Several approaches have been proposed for the
analysis of spatio-temporal processes but we prefer space-time autoregressive mov-
ing average (STARMA) models (see [7]) since these models are parsimonious and
have been widely used for the data analysis in many areas of research, such as
meteorology [9], criminology [7] and air pollution [1] etc.

The data we used in this paper is described in Sect. 3.2. Section 3.3 introduces
the multivariate regression models with STARMA errors. The model estimation,
selection of orders, and the diagnostic checking are also included in this sec-
tion. The asymptotic properties and simulations of the estimators are considered in
Sects. 3.4 and 3.5. The application of this model to the analysis of the temperatures
in the Antarctic Peninsula is considered in Sect. 3.6. The results are summarized in
Sect. 3.7.

3.2 Data

The Scientific Committee on Antarctic Research (SCAR), an inter-disciplinary com-
mittee of the International Council for Science (ICSU), established a Reference
Antarctic Data for Environmental Research project which produced a database
called READER. The database is posted on the following website: http://www.
antarctica.ac.uk/met/READER/

The database consists of monthly mean surface temperatures of several Antarc-
tic meteorological stations for recent decades. Temperatures are recorded in Celsius
scale. The monthly mean data are computed using the mean of the 6-hourly syn-
optic observations. The longitude and latitude of each station are also provided by


http://www.antarctica.ac.uk/met/READER/
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READER. The details of the database and the quality control of the observations is
discussed by [12].

There are some missing values in the database. Each missing value is estimated
by taking the average of previous two observations and next two observations in the
same month of this location. For example, the estimate of the missing value in June
1995 can be obtained by taking the average of the observations in June 1993, June
1994, June 1996 and June 1997. To obtain reliable data, we only consider continuous
periods without many missing values.

The database includes records from research stations and automatic weather sta-
tions. Since most automatic weather stations only have very short records (since
1990), we will not include these stations in our analysis. Table 3.1 shows the
location and available records of the five meteorological stations in the Antarctic
Peninsula. In our spatio-temporal analysis, we use the same period for the temper-
atures analysis at different locations. To keep the balance between longer records
and more locations, we decide to use the temperatures at Bellingshausen, Maram-
bio, Faraday/Vernadsky, Orcadas and Rothera over the period from January 1978 to
December 1998 in our analysis (the observations from January 1999 to December
2000 will be used for the prediction purpose).

Suppose we have two locations A and B, with latitude (6;, 6,) and longitude
(¢1, P2) respectively, then the great circle distance between A and B is given by:

3693.0 arccos[sin 6} sin 6, + cos ¢ cos ¢, cos(¢p; — P2)], (3.1

where 3963.0 is the radius (in miles) of the spherical earth. Therefore, the pairwise
distances of the above five stations are given in Table 3.2.

Table 3.1 The list of selected locations

No. Meteorological station Period Latitude Longitude
1 Bellingshausen Jan 1969-Dec 2008 62.2S 58.9W
2 Marambio Jan 1971-Dec 2003 64.2S 56.7TW
3 Faraday/Vernadsky Jan 1951-Dec 2006 65.4S 64.4W
4 Orcadas Jan 1904-Dec 2000 60.7S 44.TW
5 Rothera Jan 1978-Dec 2008 67.5S 68.0W

Table 3.2 Distances (in miles) between different sites at the Antarctic Peninsula

Site Bellingshausen =~ Marambio  Faraday/Vernadsky = Orcadas  Rothera
Bellingshausen 0.000 155.6 281.3 498.9 459.8
Marambio 155.6 0.000 248.3 465.5 402.1
Faraday/Vernadsky 281.3 248.3 0.000 713.7 179.3
Orcadas 498.9 465.5 713.7 0.000 860.2

Rothera 459.8 402.1 179.3 860.2 0.000
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3.3 Multivariate Regression Models with Space-Time
ARMA Errors

3.3.1 The Model

Let Y;; denote the observation at time ¢ at location i where i = 1,...,N. Let
X, = (Xi,....X,;) be a deterministic vector of covariates at time point #
t=12,....T)LetY, = (Y1s,...,Yn:) then Y, follows multivariate regres-
sion models with space-time ARMA (py,,...1,4m;...m,) €rrors if it can be written
in the form:

Y, = f(izvé) +Z,

(3.2)

P Ak O q mg o
Z, =3 2 uWOIZ, o+ 3 > 0uWWe  +g
k=11=0 k=11=0

wheret =h+1,...,T and h = max(p, q).

Though the methodology to be described here can cope with nonlinear regres-
sion model, here we will restrict ourselves to the linear regression models. For
convenience, we assume:

Xz = BX; ‘I'Zz

and we consider STARMA (pa,....4,>qmy,...m,) models for Z,.

In model (3.2), p is the autoregressive order and ¢ is the moving average order.
Ak is the spatial order of the kth autoregressive term and my is the spatial order of
the kth moving average term. The scalars ¢; and 60; are the autoregressive and
moving average parameters at temporal lag k and spatial lag I respectively. W@
is a non-zero N x N known weighting matrix at spatial lag /. Furthermore, {g,} is
assumed to be i.i.d. random vectors, with:

E(Qz) = Q
and

e, 5 =0

B = {0 20

It is further assumed that the zeros of
P Ak P Ak

det | In+ Y Y WK | and det| Iy +> > 0 wOF

k=11=0 k=11=0
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are outside the unit circle, which ensure the invertibility and the existence of a
weakly stationary solution to the space-time ARMA models given by (3.2). We also
assume that the above polynomials have no common zeros.

We have to define the weighting matrix based on the knowledge of the physical
features of the spatial locations. Since the system under study is irregularly spaced,
one reasonable approach to define the weighting matrices is to assume the spatial
orders Ay = 1 and my = 1 for all k and we have:

WO =1y and W =w

and the (i, j)th element of W is given by:

1/ l'ji?éj
» d 7 33
Wi 0 i=j (3.3)

where d;; is the distance between the sites i and j. The weights are also scaled such

N
that " w;; = 1,foreachi =1,2,...,N.
=1

Such a definition of the weighting matrix is based on a reasonable idea: the tem-
peratures from site i and j (i, j = 1,..., N) which are close to each other will be
more correlated. Hence, a greater weight of the effect of temperatures at site j will
be considered when we want to estimate the temperatures at site i.

By considering the particular weighting matrix described above and by restricting
the function f(X,, ) to be linear, the model we consider, as a special case of the

model (3.2), is given by:

Y, =BX, + 2,

p q (3.4)
Z, =) (roIn + W) Z,_ + > (Oodn +01W)e,_; + &,
k=1 =1
wheret = h+1,...,T and h = max(p,q). B is a N x r matrix of unknown
regression parameters. The By, , (im = 1,...,N;n = 1,...,r) element of the

parameter matrix B represent the effect of the nth covariate on the mth location. The
scalars ¢rg, ¢r1 and Oxg, Ok are the autoregressive and moving average parameters
at temporal lag k and spatial lag 0 and 1. W is a non-zero N x N known weighting
matrix which is defined in (3.3). {g,} is assumed to satisfy the conditions stated
earlier. To ensure that there exists a weakly stationary solution and the invertibility
is satisfied, similar conditions as model (3.2) are imposed.

3.3.2 Model Estimation and Model Selection

The model selection is done in two stages. Firstly, we choose the appropriate covari-
ates in (3.4) under the assumption that {Z,} are i.i.d. Then the residuals {Z,}
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are obtained by using least square estimation. We assume {Z .+ follows a pure
STARMA models and the autoregressive order p and the moving average order
q are determined by minimizing BIC criterion given by:

Tn|%,| 4 2k In(T) (3.5)

where k is the number of parameters in the STARMA model. T is the number of
observations and X, is the estimate of the covariance matrix of {&,} obtained by the
methods of model estimation described later.

Suppose the autoregressive order p and the moving average order ¢ have been
identified, to estimate the regression parameters By, »,(m=1,... ., N;n=1,...,r),
the autoregressive parameters ¢ro, ¢r1 (kK = 1,..., p) and the moving aver-
age parameters 09, 61 (I = 1,...,q) in model (3.4), the weighted least square
estimation procedure is used. The weighted sum of squares is defined by:

T
S= Y X lg t=h+1...T (3.6)
t=h+1

where

p q
&=Y,~BX,— > ($roIN + p1 W)X, — BX, )= > (BroIn + 01 W)e,_
k=1 =1

and X is the current estimate of the covariance matrix of &

The weighted sum of squares can be minimized by using Newton—Raphson tech-
nique. This technique provides iterative estimates of the parameters (see [6]) but
good initial estimates are still necessary. The initial estimate of regression param-
eter B is obtained by assuming that Z, is a sequence of independent random
variables. For the regression model with STAR errors, the initial estimates of autore-
gressive parameters are obtained by regressing Z, on Z,_l, WZ,_I, e ,Zt_p,
WZ t—p- For the regression model with STARMA errors, we first generate {&:}
through a regression model with high order STAR errors, then the initial esti-
mates of autoregressive and moving average parameters are given by regressing Z :
onZ, ,WZ,_,.8,_,,W_,.... The details of the procedure to find the initial
estimates for STARMA models was described in [10].

When the fitted model is obtained, it is important to test whether the residuals {&, }
are independent. The sample space-time autocovariance function of ¢, is defined by:

N

&)y
Y1k (s) = trace ([W W 28(5))

where
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The sample space-time autocorrelation function is given by:

Vik(s)

PS) = o O O

Pfeifer and Deutsch [7] suggested that specific patterns can be observed by plotting
Pro(s),(s =1,2,...) for! =0, 1, if residuals {, } are autocorrelated.

3.4 Asymptotic Properties and Simulations

Now we consider the model (3.4) described above. In this model, suppose g, =
(¢1z5-..,eNt)" and covariate vector X, = (Xi,...,Xr)'. Let w;; denote the
(i, j)th element of the weighting matrix W. The weighted sum of squares in (3.6)
is denoted by S. To derive the asymptotic properties of the least square estimates of
the model (3.4), we make the following assumptions:

e {g,} are i.i.d. with mean zero and variance 02 y.
e The zeros of

p p
det (IN + Z((bko + ¢r1 W)gk) and det (IN + Z(@ko + 61 W)gk)
k=1 k=1

are outside the unit circle.
o E(Z, 4Z, 1) isfinitefork, k' =1,..., p.

e The covariates X;; are bounded, and further for fixed i, j = 1,...,r and k,l =
1,....p
T
lim —
Tglloo Z Xi—k)X je—1)
t=h+1

exists and the r x r matrix with (i, j)th element given by lim % > XX,
T—>00 " y—pt1
is positive definite.

Furthermore, we assume X, = o021y for simplicity. Under this assumption, the
sum of squares is given by:

T
1
S=— Z gg, t=h+1...T
t=h+

Theorem 3.1. Suppose ¥ = {Bmn, Pxo, Pr1, 610,011}, withm = 1,...,N;n =
rik=1,...,p;1 =1,...,q. Theleast square estimates ¥ are asymptotically
normally distributed with mean ¥ and with covariance matrix given by:
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Cu0 0 0 o |

0 sz C23 C24 C25

0 C3; C33 C34 Css

0 Cup Cy3 Cys Cys

0 Cs; Cs3 Cs4Css

where

1 928 iy 3811
Cip= lim ———— =
N e T 0By 9B 02 P T t ;1 12; 0Bm.n 0B

withm,m' =1,...,N;n,n’ =1,...,r,and

Cor = E 2 ) —1XT:2E(Z’Z)
27 U\T dgrodgnng ) — To? G THETREH

T

1 9% 1
Cys=E[= _ BT Wz
? (T8¢k13¢k/1) To? t=2h-:|-1 (WZ,_ ) WZ, )

Crom Copm E( LS Y _ ! XT:ZE((WZ YZ, )
23 — L32 = T a¢k08¢k/1 - TUZ et Lr—k) Z—k/

withk, k' =1,..., p, and

1 %8 1 < . .
Cas=E(Fopa—]) =gz 2 2Lty +eiype)
109710 t=h+1
1 %S 1 < ) )
C55 =F 739 50, = To2 Z ZE((WQI—I) Wgt—l’ + (Wgt—l—l’) W§t)
noewr t=h+1
1 9%s ’
Cys=Css=E 73910391/1 T02 Z 2E((We,_ l) &+ We)e,)

t=h+1
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with[,l’ =1,...,q, and

1 %S [
Cyuy=Cp=E|= = 2E(Z!
= Co = E (T ) = 7o 2 M
1 s 1 <
Cis=Cs3 = E (= = S 2E(WZ, )W
35 53 (T3¢k13911) T02t=h+1 (WZ, ) Wery)

Cos=Coym E(L_OS Y ! XT: 2E(WZ,_)e,,)
25 — L52 — T a¢k03911 - TUZ et Lr—k) &r—1

CouzCporp(L 05 Y ! XT:ZE((Z Y We, ;)
TR TN T 0k 0010 T To? L TUUSTI AT

35

Proof. To prove Theorem 3.1, we follow the method of [8] given for univariate
regression models with ARMA errors. We will first show that the first order deriva-
tives of S with respect to ¥ evaluated at true values asymptotically converge to zero
and the second and third derivatives evaluated at true values converge to some con-
stants in probability. Then we verify that the estimates ¥ are consistent and evaluate

the covariance matrix by a first order Taylor expansion of the equation:

198
— =0
T gy

Finally we will establish the normality of the estimates.
As the first step to the proof for Theorem 3.1, we will show that:

—— — 0 inprobabilityas 7 — oo

where ¥ = {Bp n, bro, Px1, 010,01}, m =1,... . Nin=1,....r;k =1,...

[=1,...,q and h = max(p, q).
Differentiating S with respect of B, ,, we have:

T N

N 2 88”
=k Y Y,
0Bm.n i erwrirt 0B n

» D5
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where
»
96 Xt + Y (Prawii + dro) Xng—ky ifi=m
gir  _ k=1
0Bmn | & e
’ Y (Pawii + dro) Xng—k) ifi#m

k=1

¢j; are independent with mean zero and 3?;" L are finite and independent of ¢g,.
m.n

Consequently, from the weak law of large numbers, we have:

198
T 9B n

— 0 in probabilityas T — oo.

Differentiating S with respect of ¢y, we have:

T N

LAY 2 88”
Y Y,
ko 0% 57 15 9k
where
ae; 4
a¢: =~Yie—t)+ ) BinXna—t) = —Zig—k). forall i=1,.N.
0

n=1

N
Let M; = 0—22 > Zi—k)é&ir, using the Markov’s inequality, we have:
i=1

1 & 1 d ’
P T 2: M;| >« 58—2(T)_2E 2: M;| ., forall &> 0.
t=h+1 t=h+1

Itis easy to prove that Z; ;_x)€;; have zero mean and uncorrelated over time . Since
M; is sum of Z;_ (i)eiz, E(M;) = 0 and E(M; M) = 0 for ¢t # s and we have:

2
T T T
E{ Y M| = > EM)+2) EMM)= ) EM),
t=h+1 t=h+1 t>t’ t=h+1

for t,t'=h+1,...,T.

Furthermore, since E(M;)? < oo, we have:

|

T
zs) < SO Y EMY = ST max(EM)).

1 T
T 2 M
t=h+1 t=h-+1
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Therefore:
T
dim P | > M|ze|=0 forall £>0

t=h+1
and hence:
1 & 1 9S . .
— Z M, = — — 0 inprobabilityas T — oo, for k=1,...,p.
Tt=h+1 Ta¢k0

Differentiating S with respect of ¢, we have:

_ 381t
8¢k1 - 02 Z Z

t=h+1i=1
where

0¢eir

a¢k1 = —ZW,M(YM — Z B, an(t k)) = Zwlu u(t—k)-

u=1

Since W is a known matrix, the asymptotic properties of - 08

T 36T is similar to L 05—
Therefore, the proof will be omitted.

1
T 3¢xo
Differentiating S with respect of 6;9, we have:

0g; de;
8910 - 02 Z Z it gy where Wj; = —¢ia)

t=h+1 l—l

Since {&} is a sequence of zero mean ii.d. random variables, we have
E(gi¢-neir) =0forl =1,...,q.

We can also prove that E(g;—1&ir, &i(s—1)8is) = 0 for t # s. Following a
similar procedure described above, we can show that

——— — 0 inprobabilityas T — oo forall [ =1,...,q.
T 3659

Similarly, since we have:

0eis
89; - 02 Z Z %, eir Wwhere 89; = - Z WiuEu(t—1)-
1 t=ht1i=1 11 1

u=1
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Wwe can prove:

1 a8
——— — 0 inprobabilityas 7 — oo forall [ =1,...,q.
T 30;;

Therefore, the first derivatives of § with respect to all the parameters By, », ko,
dk1, 010, 011 converge to zero in probability.
Now we show:

1 9%s
T 0woy’

— C inprobabilityas 7T — oo

where ¥ = {Bm.n, Pko, Pk1,610. 01}, m =1,....N;n=1,....,r;k =1,..., p;
I =1,...,q and all the elements in the matrix have been defined earlier.
Obviously, we have:

1 RN

?m —> Cll in probablhty as T — oo.

For Ve > 0, by Markov’s inequality, we have:

p (| 1 RERY Cos > ) - IE (| 1 RERY C |)
S - P - e S .
T 0rodrro 2 T e T 0¢pro0¢rro 2

Since

1 RERY
T 0000

>8)=0.

C,, isdefinedas FE ( ) , we have:

1 2
lim P (‘—B—S — C22
T—o0 T d¢roddrro

Therefore, we have:

1 9°S

————— — Cy, inprobabilityas 7 — oo.
T 0¢rod¢ro

Similarly, we can prove that the other terms converge to their corresponding expec-
tations.

Differentiating S with respect to regression parameters By, , and autoregressive
parameters ¢, we have:

32—5 22 XT: ﬁ:( T + 0eir 38,’;)
8Bm,nad’ko o - 0By, na¢k0 aBm,n 8‘kaO

h+1i=1
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where

8281‘[ 88it
—— = X, 4—k) and
aBm,n 8¢k0 n= a(ka

= Zi(t—k)-

. 326‘” 381'{ : LV .
Since both B i1 and B are finite, it is easy to show that:

1 9%S

——————— — (0 inprobabilityas T — oc.
T 8Bm,n 8¢k0 P Y

Now we consider:

?s 2 XT:
aBm,na(]bkl o2 -

i ( Pei Ve asl-,)
—\0Bundprs ' B I

h+1i=1
where
828“ agil al
——— = w; X,,¢u—k) and = WinZy(t—k
0B11,0 0¢r1 n= 01 ; =0
and w;; is the (i, j)th element of weighting matrix W for (i,j = 1,...,m).
Obviously, the asymptotic properties for 33;32% is similar to zm,?,z,,—sétme» SO we
have:
1 928
————— — 0 inprobabilityas 7 — oo.
T 3Bm,n 0Px1
Since:
928 2 & 0%ei; dej; 0Ogis
R A0 2 Z Z it + e
0B 000 o ‘ 0B, 06010 0B 39;0
’ t=h+1i=1 ’ ,
where
8281‘[ 881‘[
——— =0 and — = —¢g;y—
9Bm.n 010 30 He=h

by using the weak law of large numbers, we can easily show:

1 928 1 9S

=7 —0 d =———F+——0 i babilit T .
T 3B 310 — an T 3B 900 — 0 in probability as — 00
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Therefore, all the second order derivatives converges to matrix C which is given
earlier.

Similarly, it is not difficult to prove that the third order derivatives of S are
uniformly bounded, i.e., we have:

! ) M i babilit
——————— — M, i, inprobabili
T 0w, 0w, 0w, o oves 1P Y
where ¥ = {Bmn, dr0,Pr1,00,.61}, m = 1,...,N;n = 1,...,r;k =
IL...,p;l=1,...,qand iy, iz, iz =1,...,mn + 2(p + q). M;,i,i; is a constant
independent of 7.

Now we derive the covariance matrix of the estimators.
_ For convenience, let u =mn + 2(p + ¢). Suppose ¥ is the true parameter and
¥ is the least square estimate, also ¥* lie close to the true parameter. The Taylor
expansion for the first order derivatives of S with respect to ¥ is:

T35 b= 775 +( A B LY A
Tow V=" T g YTV owaw Y= ToT ' W(?;)
where the i;th row of Q is given by:
RERY
U -y, - -
( 0) T 0W;, 09,09,

We have shown earlier that the first order derivatives on the right hand side of
(3.7) converge to zero in probability and the second and third order derivatives con-
verge to constants. Hence, by a theorem of [3], we have a consistent least square
estimate of ¥ with probability approaching 1 as T — co. Because the consistency
of ¥, 0 in (3.7) converges to zero. We also know that:

1aS 1 3*S
T3w lg—g=10 and T 3030 |g—g,— C in probabilityas 7T — oc.

Hence, from (3.7), we have:

1 aS R
ﬁﬁ lo=w, +«/T(l1/ —Y)C—>0 as T — oo.
Therefore:
1,08 N C e
ﬁC P2 lg—w, and T (¥ —Y¥%) have the same asymptotic distributions.

Since we have shown earlier that the expectation of % g—i |w=w, equals zero, (lf/— )
will also have asymptotically zero mean. The sample covariance matrix of ¥ is
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given by:

_ 10SaSY\ .,
=—=C im -
T T—o00 T 0¥ 0¥

It is straightforward to show that:

105 9dS 1 028
E (Tﬁﬁ) =25 spaer)

this implies:
1 2
r=_chwc!=_ch
T T

We now establish the asymptotic normality of the estimates. We will first study
the asymptotic normality of the regression estimates B,,,(m = 1,...,N;n =
1,...,r). We note that ai’;’ﬂ are finite and independent of ¢&,. Therefore, 3 gS
is the sum of independent random variables with different variances. Since we have
, it follows that:

ﬁmte

881'[ 881[
1r5nza§XT (3Bm,n) Z Z( ) —0 as T — oo.

t=h+1i=1

Hence, the Lindeberg condition is satisfied and \/LTaBa% are asymptotically nor-

mal by using the Lindeberg Feller central limit theorem. Since J;TI -1 3 gms.n and

VT (B — Bgn.nyo) (Where Bpnyo is the true value of the regression parameter)
have the same asymptotic distribution, it is straightforward to conclude that the esti-
mate of regression parameters By, 5 is also asymptotically normally distributed and
any linear combination % > em naBa—S is also normally distributed. Therefore,

\/L a5 1s distributed asymptotically as a multivariate normal.

We now establish the asymptotic normality of the autoregressive and moving

average estimates. From the previous proof, all the terms a‘;‘z 5 32&1 , 33050 880% are

linear sums of stationary and uncorrelated series. Moreover, they are linear sums of
: : 1 1 a8 1 9S8
martingale differences. Hence, by a theorem of Billingsley [2], T 360" JT

\IF 33050 ‘/L 36, are asymptotically normal. Since any linear combination of above

terms are asymptotically normal, the first order derivatives of S with respect to
the autoregressive and moving average parameters are asymptotically multivariate
normal.

Therefore, the least square estimators of all the parameters are asymptotically
multivariate normally distributed and hence the results of Theorem 3.1. O
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3.5 Simulation Study

The estimation methodology described above is used for simulated data. All the sim-
ulations were performed using pseudo-normal random numbers. Space-time data
sets were simulated from different regression models with different STAR(1;) and
STARMA(1y, 1) errors. We consider a system of five locations with the weighting
matrix given by:

0.0000
0.4260
0.2441
0.2984
0.1907

0.4539
0.0000
0.2766
0.3199
0.2181

0.2510
0.2669
0.0000
0.2086
0.4892

0.1415
0.1424
0.0962
0.0000
0.1019

0.1536
0.1648
0.3831
0.1731
0.0000

This weighting matrix is calculated based on the real locations of the five sta-
tions (Bellingshausen, Marambio, Faraday/Vernadsky, Orcadas and Rothera) in the
Antarctic Peninsula.

3.5.1 Model 1

We have generated space-time data set from the model given by (the number of
locations is assumed to be 5):

Y, =BX, + 2,

~ MN(0, I
Z,=(04Iy —02W)Z,_; +¢, & (0.15)

(3.8)

where:
3.9)

and

2.0
1.5
2.0
1.5
2.0

3.0
3.0
3.0
3.0
3.0

3.0
3.0
3.0
3.0
3.0

B=| -50
5.0

5.0

(3.10)

3.5.2 Model 2

We have also generated another time series from a regression model with STARMA
errors which is given below:
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Y, =BX, + 2,
Z,=(04Iy -02W)Z,_, t=2,....T & ~MNQ,Is) (3.11)
+(03Iy +02W)e,_ + &

where the covariates and the regression parameters B are same as in model 1.

3.5.3 Model 3

Besides, we have generated time series from the following regression model with
STARMA errors:

Y, =BX, + 2,
Z,= 03Iy +01W)Z,_, t=2,....,T & ~MNQ,Is) (3.12)
+(02Iy —01W)e,_; + ¢,

where:

t 2xt . 2wt
X, =11, =, cos —,sin —
T 12 12

and

=50 2.5 3.0 30
=50 2.5 3.0 30
B=] -50 25 30 3.0
=50 2.5 3.0 3.0
=50 2.5 3.0 30

Tables 3.3, 3.4 and 3.5 show the average estimates of the parameters of interest
from 100 replications for each T in the above three models. From the results of
the tables we observe that the accuracy of the estimates increase with the sample
size for all the models. Moreover, the standard deviations (given in the bracket of
estimates) of all the estimates, as expected, decrease with the sample size. Therefore,
the simulation results shows that the estimates are asymptotically consistent.

3.6 Real Data Analysis

In this section, we consider the analysis of the temperatures of Antarctic Penin-
sula by using multivariate regression models with space-time ARMA errors. The
data described earlier consists of 252 monthly mean surface temperatures recorded
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Table 3.3 Simulation results for model 1

X. Gao and T.S. Rao

Sample size T =100 T =200 T = 400
B 2 1.9998(0.1605) 2.0064(0.1224) 2.0047(0.0872)
]§2_2 1.5067(0.1948) 1.4951(0.1157) 1.5061(0.0934)
]§3_2 2.0317(0.1917) 1.9782(0.1200) 2.0039(0.0859)
]§4_2 1.4873(0.1724) 1.4825(0.1170) 1.4977(0.0854)
]§5_2 1.9980(0.1795) 2.0137(0.1150) 2.0122(0.0947)
qfvm 0.3969(0.0114) 0.3978(0.0084) 0.3994(0.0055)
q311 —0.2021(0.0207) —0.1991(0.0150) —0.2022(0.0113)
Table 3.4 Simulation results for model 2

Sample size T =100 T =200 T = 400
B 2 2.0159(0.2168) 2.0067(0.1472) 2.0044(0.1022)
éz.z 1.5161(0.2238) 1.5122(0.1801) 1.4990(0.1086)
é}_z 2.0042(0.2458) 2.0032(0.1552) 2.0067(0.1099)
I§4_2 1.5133(0.2016) 1.5013(0.1535) 1.4910(0.1005)
]§5_2 2.0233(0.2467) 1.9952(0.1604) 1.9822(0.1102)
qAﬁlo 0.3963(0.0203) 0.3966(0.0144) 0.3974(0.0095)
q31 1 —0.2027(0.0370) —0.2020(0.0293) —0.1997(0.0183)
ém 0.3016(0.0223) 0.3028(0.0148) 0.3024(0.0091)
él | 0.2035(0.0363) 0.2014(0.0255) 0.2005(0.0157)
Table 3.5 Simulation results for model 3

Sample size T =100 T =200 T = 400
B 2 2.4904(0.1832) 2.4979(0.1093) 2.4938(0.0723)
éz_z 2.5080(0.1694) 2.5050(0.1363) 2.4947(0.0792)
é}_z 2.5066(0.1665) 2.4788(0.1240) 2.4965(0.0842)
I§4.2 2.5143(0.1731) 2.4915(0.1187) 2.5049(0.0792)
]§5_2 2.5015(0.1677) 2.5106(0.1440) 2.4969(0.0811)
qAﬁlo 0.2961(0.0268) 0.2989(0.0207 ) 0.2974(0.0146)
q311 0.0995(0.0451) 0.0997(0.0330) 0.1062(0.0228)
ém 0.1990(0.0274) 0.1990(0.0209) 0.2020(0.0159)
él | —0.0994(0.0489) —0.0983(0.0365) —0.1043(0.0253)

from January 1978 to December 1998 at the five meteorological stations (Belling-
shausen, Marambio, Faraday/Vernadsky, Orcadas and Rothera) The time series plots
of the temperatures for each station is given in Fig. 3.1. All the plots suggest strong
seasonality of the series with period 12 months.

It is reasonable to use the inverse distance between two sites as the correspond-
ing element of the weighting matrix since the correlation between the residuals

{Z,} after fitting the linear regression model in (3.13) from two sites seems to be
inversely related to the distances (see Fig. 3.2). The corresponding stations of each
correlation are shown in the label of each point and the number of each station
is given in Table 3.1. For example, the point labeled (2, 4) represents the distance
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Fig. 3.2 Correlation between two series and the corresponding distance of the two sites
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between Marambio and Orcadas and also the correlation between the residuals from
the above two sites after fitting linear regression models.

The weighting matrix is calculated by the Definition 3.3, then it is standardized so
that all rows add up to one, thus the weighting matrix W given in (3.8) is obtained.
We use the following multivariate regression model with space-time ARMA errors
considered earlier to fit the data at the five locations of Antarctic Peninsula:

Xt = BX; +Zt

P q 3.13
Z, = 3 Groly + $aW)Zyp + 3 Groly + 0nW)e, , +e, 1
k=1 =1

where Bsx4 is a matrix of parameters that need to be estimated. We have:
E (Qz) =0

and

E(giglys) = 02’8’ s £ 8

Since the temperature series of all the five locations have mean and trend and
moreover they show strong seasonality with period 12, we use covariate vector
X, =11, %, cos %, sin % ’. Other assumptions of this model are same as (3.4).
The autoregressive order p and the moving average order g are determined by min-
imizing BIC criterion. From the results in Table 3.6, we observe that the BIC is
minimum when p = land g = 1.

By using the estimation procedure described before, the estimates and corre-
sponding standard errors (shown in the bracket) are calculated and are given in the
following matrix:

—2.4882(0.1723) 0.3418(0.2925) 3.1384(0.0829) 2.6238(0.0835)
—8.5167(0.2403) 0.0646(0.4089) 5.6156(0.1267) 2.2886(0.1276)
B = | —4.5733(0.1807) 2.2394(0.3070) 3.0881(0.0896) 3.5517(0.0900)
—3.6615(0.1875) 0.5901(0.3186) 4.2916(0.0943) 3.1598(0.0949)
—5.7757(0.1978) 2.2595(0.3362) 4.4711(0.0993) 4.0392(0.3169)

Table 3.6 Model selection for STARMA models

Order p=20 1 2 3

q=0 705.9 715.8 724.2
1 695.7 667.2 678.2 684.5
2 679.5 674.9 680.7 681.3

3 686.5 685.2 680.7 691.3
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Fig. 3.3 Space time ACF for residuals {2, }

$10 = 0.3374(0.0607) 1 = 0.2594(0.0727)
f10 = —0.0119(0.0541) 617 = —0.1084(0.0559).

The results in the above table show that the estimates of mean (first column
of E) and harmonic components (third and fourth column of B) at all the loca-
tions are significant. The trends (second column of B) are significant at level 1%
at Faraday/Vernadsky and Rothera. The trends are also significant at level 10% at
Orcadas but they are not significant at other locations. This analysis confirms that
the warming at Faraday/Vernadsky and Rothera are very significant, compared to
other stations. The results also show that the estimates of autoregressive parameters
and the moving average parameter at spatial lag 1 (éll) are significant.

Figure 3.3 shows that the residuals {é;} obtained from the fitted model are
not autocorrelated since autocorrelations at most lags are not significant and there
are reasons that they are normal and hence the assumption that the residuals are
independent is satisfied.

After fitting the model to our temperature data, we are interested in forecasting
the temperatures of each site in the future. Let Y, be an vector of observations at
each site at time . Let Y, (/) be the predictor of Y, ; made at time ¢. The mean
square error of the / step predictor is given by:
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-12
0
Fig. 3.4 Predictions of the temperatures from January 1999 to December 2000

M) = E[X.()-Y, )]

The optimum predictor of Y, , ; which minimizes the mean square error is:

~

L-‘rl = E(ZI+Z|L’LL‘—19"~-)~

For our model, the one step predictor for the temperatures is given by:

Yii1 = BXi1 + ($rols + puuW) (Y, — BX,) + (Brols — 911W)§t-
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The one step prediction for the temperatures from January 1999 to December
2000 at the five stations are compared with the real data over the same period. The
comparisons of the predictions with the true values are given in Fig. 3.4 (the dash
line is the prediction line). The results show that all the predictions are close to the
true observations.

3.7 Conclusion

In this paper we examined the temporal and spatial variability of the temperatures
at the five locations in the Antarctic Peninsula. Besides fitting linear regression
models to temperatures, we consider multivariate regression models with space-
time ARMA errors to study the spatio-temporal variation. From our analysis, the
monthly mean surface temperatures at all the locations in the Antarctic Peninsula
are increasing over the period from January 1978 to December 1998. The largest
warming trends are found at the Faraday/Vernadsky and Rothera, with the warming
rate of 1.0664 °C/decade and 1.0759 °C/decade respectively. The trends at the above
two locations are also significant at 1% level. The temperatures at Orcadas is also
increasing at 10% significance level, with the warming rate of 0.5901 °C/decade.

A warming rate of 1.01 °C/decade at Rothera over the period from 1978 to 2000
is also reported by [12]. However, the estimated trend given in [12] is not signifi-
cant because of the lack of efficiency by using simple linear regression models to fit
temperature data. From our analysis, we believe the substantial warming occurred
at Rothera station is significant at level 1% since the seasonality and temporal
autocorrelations have been taken into account.
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Chapter 4
The Generalized von Mises—Fisher Distribution

Riccardo Gatto

Abstract In this chapter we introduce the broad class of generalized von Mises—
Fisher (GVMF) distributions on the unit hypersphere S”~!, which arises from a
generalization of the von Mises—Fisher distribution. This class of distributions has
some important information theoretic properties. It is shown that, under constraints
on some moments along some fixed directions, and using Kullback—Leibler infor-
mation as measure, the closest distribution to any predetermined one on S?~!, has
the GVMF form. Lower bounds for the Kullback-Leibler information in this con-
text are also provided. Several connections between GvMF and other directional or
linear distributions are given. GvMF distributions can be re-expressed in terms of
generalized von Mises distributions when p = 2. GvMF distributions arise as offset
multivariate normal distributions, as offset singular normal distributions and as off-
set distributions from an exponential spherical type of distributions. Various forms
of GvMF densities which feature uni- and multimodality, with different shape of
modes, girdle and with other particularities are graphically illustrated.

4.1 Introduction

In this chapter we introduce the generalized von Mises—Fisher (GvMF) direc-
tional probability distribution, motivated from its high flexibility, its information
theoretic properties and its characterizations in terms of other important distri-
butions. Directional data which are directions in R?, p = 2,3,..., and can be
represented by points on the surface of the (p — 1)-dimensional unit hypersphere
SP~1 = {x € RP|x"x = 1}. Most practical situations are for circular data, where
p = 2, and for spherical data, where p = 3. Many results of this chapter are given
for the general case of p > 3, because it is conceptually similar to the case of p = 3.
Spherical data arise in various fields such as physics, astronomy and earth sciences
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and can have experimental or observational origins. A probability distribution on
SP~1 is a directional probability distribution. Two general references on directional
statistics are [10] and [6]. In the following we are only concerned with absolutely
continuous directional distributions with respect to (w.r.t.) the uniform distribution
on S?~1. When p = 2 it is generally convenient to represent a random unit vector
by its angle w.r.t. a chosen null direction and with the radian measure, for example.
This random angle has a circular density, w.r.t. the Lebesgue measure on S, which
is a non-negative 2x-periodic function on R integrating to one on [0, 27). In gen-
eral we can re-express x € R?\{0} in angular coordinates by the diffeomorphism
x = g(r,01,...,0,-1) which, for p > 3, is given by

x1 =rsinfysinb,...sinf,_»cosf,_1,

Xy =rsinfysinb,...sin6,_5sinb,_q,

Xp—1 = rsinf; sin by,

Xp = rcosby,

wherer > 0,0 < 60; <mfori =1,...,p—2and0 < 0,_; <2n.Forp = 3,0, is
the co-latitude and 65 is the longitude. The determinant of the Jacobian of g is given
by J(r;61,...,0p—2) = Pl ]_[f;; sin?~/ 0;_1,for p > 3, and it is equal to r for
p = 2. Jacobi’s theorem yields that for any continuous function f : SP~! — R,

[, fedow -
2m T 2w
/ / f(g(l;(?l,...,(9,,_1))J(1;(91,...,9p_2)d(91...d@I,_I,
0 0 0

where do (x) denotes the infinitesimal surface area on S?~! and around x on S?~1.
From this, the surface area of S?~!isa, = Jsp—1do(x) = 2P/ (p/2), T
denoting the gamma function. Hence, the uniform or isotropic probability distri-
bution over P~ is U[A] = [, do(x)/ap, for A C SP~1 which is the unique
invariant distribution under rotation and reflection. For a probability distribution P
over S”~! and for A C SP7!, P[A] = [,(dP)/(dU)dU, where (dP)/(dU) is
the Radon—-Nikodym derivative or density of P w.r.t. U. All directional densities
presented in this chapter are given w.r.t. U.

We now introduce a class of probability distributions on S?~! which generalizes
and unifies several other existing distributions. The generalized von Mises-Fisher
distribution of order k (GvMFy,) is the probability distribution P on S?~! having
the density w.r.t. the isotropic distribution U at x € S?~!

def dP

xX|lmq,....my;c1, ... Chilit,...,if) = —
Sf(x|m, my; ¢y ki1 k) J0

k
(x) o< exp g > Cr(miz)"’§ :

r=1

“.1)
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where m, € SP71, ¢, e R, forr = 1,...,k, iy < ... <ix € {1,2,...} and, to
avoid over indentification, 1, (mx)'!, ..., (m; x)'* are linearly independent func-
tions of x € S?~!. We denote as GVMF,(my,....my;c1,...,Cksit,...,0k) a

representative random direction of R? with density (4.1). The particular case of
k = 1andi; = 1in (4.1) yields the von Mises—Fisher (vMF) density which plays
an important role in directional statistics (see, e.g., [10, p. 168]), and we denote as
VMF(m,;c1), m; € R?, ¢y > 0, a representative random direction of R? with this
density. Thus, a GvMFy with iy, ..., known belongs to the most general expo-
nential directional distribution of [2], with density proportional to the exponential
of polynomials in x € S?7!. The reparametrization (4.1) leads however to intu-
itive interpretations of the parameters, to information theoretic optimalities and to
various characterization properties in terms of other important distributions.

In Sect. 4.2, we show that GVvMF distributions possess some important informa-
tion theoretic properties. We first show that for some fixed moments along some
predetermined directions, the closest density, in the Kullback—Leibler sense, to a
given density fj, all of them on S?~!, has the GYMF form (4.1). We also provide a
lower bound for the Kullback—Leibler information under these moment restrictions
as well as the entropy of GvMF distributions. In Sect.4.3, we show that GVYMF
distributions are closely related with other important directional and linear distri-
butions. When p = 2, any GvMF distribution admits a generalized von Mises
(GvM) reparametrization. The GvM distribution was introduced by [5]. GvMF,
distributions are closely related to the Fisher—Watson and the Fisher—Bingham direc-
tional distributions. GvMF distributions arise also as conditional offset distributions
from the multivariate normal, the singular normal and from a type of spherical
exponential distributions. Still in Sect. 4.3, we show various graphs of GvMF densi-
ties, which feature uni- and multimodality, different modal shapes, girdle and other
interesting forms.

4.2 Information Theoretic Results for Directional
Distributions

In this section we derive some optimal directional distributions w.r.t. directional
versions of Kullback-Leibler information and entropy, respectively defined by (4.2)
and (4.3) below. Under both criteria, either GVMF distributions or other directional
distributions having a GvMF part turn out to be the optima. An important con-
sequence is that these distributions are optimal solutions of a constrained prior
selection problem of Bayesian statistics. In our setting, an optimal directional prior
distribution is obtained by maximizing the entropy given some prior knowledge on
the moments along some directions.

The Kullback-Leibler information was introduced by [8] and plays a central role
in information theory. We suggest defining this quantity on S?~! also. Suppose P
and Q are two probability measures on S?~! w.r.t. the uniform distribution U and
that P is absolutely continuous w.r.t. Q, which is denoted P < Q. Then we define
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_ a S(x)
I(P|Q) = /;p_l log EdP = [Sp_l log @f@)dU(Q, 4.2)

where f and g are the directional densities of P and Q respectively w.r.t. U and
where 0log 0 = 0 by assumption. /(P|Q) is the mean logarithmic likelihood ratio
w.r.t. P or mean information per observation of P for discriminating of Q from P.
The Gibbs inequality implies that /(P |Q) is positive semi-definite, i.e.,

1(P]Q) =0, (4.3)

for all P and Q such that P <« Q, with equality iff P = Q as. I(P|Q) is
sometimes referred to as relative entropy or Kullback-Leibler distance, even though
I(P|Q) is not a metric: it violates the symmetry and the triangle rules. A related
quantity is the entropy of [12], which we suggest extending to S?~! by

dP
1)y == [ woefgar == [ g ) f@dU)

where 0log0 = 0 is assumed. H(P) measures the uncertainty inherent in P or
in f. Equivalently, H(P) measures the expected amount of information gained on
obtaining a direction from P, based on the principle that the rarer an event, the more
informative its occurrence. We denote alternatively I( f|g) for I(P|Q) and H(f)
for H(P).

We denote by DM, the rth directional moment condition on the density f given
by

[, v = o,

wherei; <...<ip e{l,2,...},m, € SP7l g, eRandr =1,...,k.
All densities in the next theorem and corollaries are on S?~! and w.rt. the
isotropic distribution U on S7~!.

Theorem 4.1. (a) The directional density [ which satisfies DMy, forr = 1,...,k,
and which minimizes the Kullback—Leibler information 1(f|fo), w.rt. a given
directional density fy, is proportional to

k
exp { > e (mﬁ&)i’} fo), (4.4)
r=1

forx € Sp-1 The parameters cq, . . ., Ck are the solutions in vy, ..., Vg of

a0
F KWi,...,vismy, .. ,mysin, .. ik fo) =ar, r=1,...,k, 4.5)
Vr

where K = log M and
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k

M@y, .. ovesmy, . myin, kS fo)=/S lengz vr(miz)i’§fo(£)dU(£).
.

r=1
(4.6)
(b) For any directional density f satisfying DM,, forr = 1,...,k, we have the
following lower bound on the Kullback—Leibler information,

k
I(f1fo) = —K(cr,....ciimy....ompiiv....igs fo) + Y cjoy. (A7)
j=1

with equality iff f is proportional to (4.4).

Proof. The basic idea of th_is proof'is due to [7].
(a) Define t,(x) = (m,x)",r = 1,...,k. In order to minimize /(f|fo) w.r.t. f
and under DM,, forr = 1,...,k, we minimize w.r.t. f the Lagrangian

k
Ln= [ glog R —Vo} SU)

= [ v hwave.,

where

k
v(g) = (logq = vty — Vo) q

r=1

and ¢ = f/ fo. Define

k
qozexp§Zvrtr+vo—1}.

r=1

We note that ¥(q0) = —qo. ¥'(q) = logq — Yk, vetr —vo + 1, ¥/(qo) = 0,
¥"(q) = 1/q. For any function 1 on S?~! between g and go, we have

L= [ [pa0) + 19w - g0@iv o)
+ 240~ 400" () | /o) U )

[ @ h@aUw+3 [ a0 - 0w W@ AWdU.
Ssr—l1 sp—1
(4.8)
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This implies

k
Lnz= [ ew { > vrte(@) +vo - 1} fo@dU),  “9)

r=1

with equality iff the second integral in (4.8) vanishes, which happens iff

k
f= exp{Zvrtr + v — 1} fo-

r=1

From the definition (4.6) and because f integrates to one w.r.t. U on S?~!, we have
et = MY(vi, ... viimy. .o mycin, ks fo). Hence we obtain

k
fx) = M_I(Vl,---,vk;ml,---,mk;il,m,ik:fo)eXP{ZVrlr(l)} Jo(x).
r=1

(4.10)
From DM,, r = 1,...,k, follows that cy, ..., cg are the solutions in vy, ..., vg of
4.5).
(b) From (4.8) and (4.9), we obtain that for any density f satisfying DM,, r =
1,...,k, (4.7) must hold and it must hold with an equality iff we take the optimal
f given by (4.10). O

Corollary 4.1. (a) For any k € {1,2,...}, the directional density [ maximiz-
ing the entropy H(f) under DM,, r = 1,....k, is the GYMFy(m,, ..., my;

Cly... Ckii1,...,ir) density. The parameters cy,...,cr are the solutions in
Vi, ..., Vg Of
a . o q 2 .
WK(vl,...,vk,ml,...,mk,tl,...,lk,1)=otr, 4.11)
r
r=1,...,k.

(b) If f is a directional density satisfying DM, r = 1,...,k, then

k
H(f) S K(c17'-‘7ck;m]7'-‘7mk;i1""aik;1)_Zcrar’
r=1

with equality iff f is the GVWMFy(my, ... ,my;c1,...,Ck;i1, ..., i) density.

Proof. (a) By choosing fo = 1 a.s.on SP~!, we find I(f|fo) = —H(f). Hence
maximizing H(f) w.r.t. f under DM,, r = 1,...,k, is equivalent to minimizing
I(f|fo) w.rt. f under these constraints. It follows from Theorem 4.1(a) that this
minimum is attained when f is a GvMFy density. The parameters cy, . . ., ¢y Which
satisfy the constraints are given by (4.11), which is exactly (4.5) with f, = 1.

(b) This part follows directly from Theorem 4.1(b). O
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As already mentioned, Corollary 4.1 can be used for prior selection in Bayesian
statistics. An appealing property of the GvMF class of distributions is its closure
w.r.t. directional moment restrictions in the following sense. All densities are again
given w.r.t. the isotropic measure U on S?~!. The proof of the following corollary
is direct and omitted. In the next corollary, we relax the inessential restriction on the
orderingiy < ... <i; <ij41 <...<ij4k.

Corollary 4.2. (a) Denote by fo the GYMF;(my,... ,m;;c1,...,cjiit, ... i)
density. Then the closest density f to fy, in the sense of minimizing 1(f| fo),
which satisfies DMy, r = j + 1,...,j + k, is the GYMF; yx(my, ... . m; x;

Clye s Cjtkiils ..., 1j+k) density. The parameters cji1,...,Cj4+k are solutions
iNVjt1, ..., Vjyk of
WK(Vj+17"'7Vj+k;mj+1’"'amj+k;ij+l7'-~7ij+k;f0) = Uy,
-

r=j+1,...,j +k.
(b) For any density f satisfying DM,, r = j +1,...,] +k,

j+k
I(f 1) ==K (Cjgte o Cipki Mgyt O+ D Cr,
r=j+1

with equality iff f is the GYMF . density given in (a).

4.3 Other Properties

In this section we first provide some other properties of GvMF distributions, which
concern the invariance, the clustering around the mode and the antipodal symme-
try, and we show a few of the various possible shapes taken by GvMF densities.
Then we show various relationships between GvMF and other directional or linear
distributions.

The first property of GvMF distributions is their invariance under orthogo-
nal transformations. If X ~ GvMFg(m,,....my;c1,...,cksi1,...,0ix) on §p1
and A4 is a p x p orthogonal matrix, then ¥ = AX ~GvMFy(Am,,..., Amy;
Cly...,Ck;i1,...,ir), meaning that the class of GvMF distributions with fixed
C1s...,Cgand iy, ..., I, is invariant under the group of orthogonal transformations
on R?. As practical consequence, the choice of coordinate system is irrelevant.

Regarding the shapes taken by the GVMF densities we can provide the follow-
ing general statements. If i, is even, then the clustering around both directions
m, and —m, increases as ¢, increases, r = 1,...,k. If iy,...,i; are even, then
(dP)/(dU)(x) = (dP)/(dU)(—x) Vx € SP~!, meaning that we have invariance
under x —> —x,x € § =1l e antipodal symmetry, and we have thus a distri-
bution for axial data. In general, the larger the values of ¢y, ..., ¢k, the larger the
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Fig. 4.1 GvMF,((0,0, 1);
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clustering around m, ... ,m;. lf my = ... = my and cq,...,cx > 0, then the
maximum of the density is at m; and the minimum at —m ;. If m;, = ... = my,
then we have the rotationally symmetry around m,. Clearly, the GvMF distribu-
tion with ¢y = ... = ¢x = 0 is the isotropic distribution. Any GvMF;, distribution
withi; = ... = ix = 1 is simply the vMF(||v]|~v; ||v||) distribution, whenever
v=Yr_ cm, #0.

Figures 4.1-4.12 show some particular GYMF densities on S2. The densities
are plotted around the unit sphere and up to unspecified proportionality constants.
GvMF, densities with i; = 1,3 and 7 are respectively plotted in Figs. 4.1, 4.2 and
4.3, all other parameters being held fixed. These figures show hence the effect of
the increase of i1 in the GVMF; density. Figures 4.2 and 4.4 have GvMF; densities
with ¢; = 1 and 2 respectively, all other parameters being equal. Figures 4.4 and
4.5 are GvMF] densities with i; = 3 and 4 respectively, all other parameters being
equal. Both Figs. 4.5 and 4.11 show antipodal symmetries, yielding distributions for
axial data. Figure 4.5 has i; = 4 even and has hence a mode and a similar antimode.
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Fig. 4.3 GvMF,((0,0, 1); i -
157) 15 ..o o
14. ...
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Fig. 44 GvMF((0,0,1); ,
2;3) Sy

Figure 4.11 is a GvMF3 density with i; = i, = i3 = 4 and hence with three
modes and three similar antimodes. Due to the choice of the other parameters, all
six modes are in fact similar. Figures 4.6 and 4.7 show two GvMF, densities and
Figs. 4.8-4.11 show four GvMFj3 densities. Finally, Fig. 4.12 shows a girdle shaped
GvMF, density.

Another existing generalization of the GvMF distribution is the Fisher—-Bingham
distribution, whose density at x € S?~! is proportional to

exp{em'x + x"Mx},

where ¢ > 0,m € SP~! and M is a p x p symmetric matrix standardized to have
null trace (see, e.g., [10, p. 174]). The restriction to matrices with null trace affects
the normalizing constant only. If we restrict M to have rank one and relax the null
trace condition, then both symmetry and unitary rank conditions are equivalent to
the existence of a vector ¢ € R” such that M = aa’. By setting a = ,/cam,,
where ¢, > 0 and ||m,|| = 1, we see that the GVMF, distribution with i; = 1 and
ip = 2 is the special case of the Fisher—Bingham distribution with rank{M} = 1.
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Fig. 4.5 GvMF,((0, 0, 1);
2;4)

Fig. 4.6 GVMF,((0, 1,0).
(0,0,1);1,1;3,5)

This latter GvMF, distribution is in fact known as Fisher—Watson distribution (see
[10, p. 177]).

Fig. 4.7 GVMF,((0, 1,0).
0,0,1);1,1;3,10)
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Fig. 4.8 GvMF;((1,0,0).
(0,1,0),(0,0,1);1,1,1;3.3,3)
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Fig. 4.9 GVMF;((1,0,0).
0,1,0),(0,0,1); 1, 1,15 15,15, 15)

For p = 2, a broad class of circular distributions which plays an important role
w.r.t. the information theoretic quantities defined in Sect. 4.2 is the generalized von
Mises of order kK (GvMg). In terms of the angular coordinate and w.r.t. isotropic or
the Lebesgue measure on S!, the GvMy, density is given by

f(9 | HUiseeos Ri3KT, oo KE) =
1 k
exp Kkpcosr( —up)e . (4.12)
271G((,k)(81,...,Sk_l;/q,...,/ck) r;

for 6 € [0,2x), kr > 0, uy €[0,21t/r),forr = 1,...,k, and where
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Fig. 4.10 GvMF;((1,0,0),
(0,1,0), (0,0, 1); 1, 1, 1; 15, 15, 15)

Fig. 4.11 GvMF;((0,0, 1),
0,1,0),(0,0,1);2,2,2;4,4,4)

Fig. 4.12 GvMF,((0, 1,0),

(1,1,0)/4/2,(1,0,0), (1, =1,0)//2;
1,1,1,1;6,6,6,6)
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k
GG ik k) =
1 2

T expi{ky cos 6 + kpcos2(0 4+ 81) + ... + kp cosk(0 + Sx—1)}d0,
T Jo

with §; = (1 —p2)modm, 8, = (w1 —pu3)mod(27/3),...,8k—1 = (1 —ur)mod
(27/ k). We denote by GvMy (i1, ..., ik: K1, ..., K ) a Tepresentative angle with
the density (4.12). This class of distributions was introduced by [9] and [5] as a
generalization of the circular normal or von Mises (vM) distribution, whose density
is given by

FO 1) = expik cos(6 — )},

1
21y (K)
for 0, € [0,2m), k > 0, where I,(z) = (2m)~! fozn cosnb exp{zcos0}do, z €
C, is the modified Bessel function of the first kind and integer order n (see, e.g.,
[1, p. 376]). Compared to the vM, which is circular symmetric and unimodal, the
GvM; distribution allows already for higher flexibility in terms of asymmetry and
bimodality.

When p = 2, many GvMF;, densities with are indeed GvMy, densities. The
following result we illustrates this fact for the case k = 3 andi, =r,r =1,2,3.

Result 4.1. Consider X ~ GvMFz(m;,m,,m5;c1,¢2,¢3;1,2,3) on S and define
6 = arg{X}. Then 8 ~ GvM3(u1, 2, 13; k1, k2, k3), where for v, = arg{m,},
r =1,2,3, and arctan : R — (—n/2, 7/2), we have

4cq sinvy 3c38inv
u1= | arctan ! 1+ 96 3 +ml{4cy cosvy + 3c3cosvz < 0} | mod 2,
4cqcosvy + 3c3cos v
(4.13)

K1 = {c? + (9/16)c§ + (3/2)cic3 cos(vy — v3)}%, (4.14)

U2 = Va,k3 = c2/2, 03 = v3 and k3 = c3/4.

Proof. Let us define 6 = arg{x}, where x € S!, v, = arg{m,} and 6, = 6 — v,
for r = 1,2,3. From mix = cos 6y, (mhx)* = cos*6, = (1/2)cos26, + 1/2
and (m}x)*® = cos® 03 = (1/4)cos363 + (3/4) cos B3 and from the fact that the
transformation from Cartesian to polar coordinates has Jacobian one, it follows from
(4.1) that the density of arg{ X} at 6 is proportional to

3 1 1
exp {cl cos(8 —vy) + ZC3 cos(6 —v3) + 562 cos2(6—vy) + ZC3 cos 3(9—1}3)}.
(4.15)
The first two summands of the above exponent can be re-written as

3 . . 3.
cosf | cicosvy + ZC3 cosvs ) +sinf | ¢y sinvy + ZC3 sin v3

= cos 6 (k1 cos 1) + sinf (kqsinpg) = xqcos(6 — py),
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with @1 and «7 given by (4.13) and (4.14). From this follows the GvM3 density at 6

1
270G (81, 82: k1, k2, k3)

expi{xy cos(0—p1)+ro cos2(0—pa)+k3 cos3(0—us3)},

where w1 is given by (4.13), k1 is given by (4.14), uo = va,kx = ¢2/2, 43 =
v3,k3 = ¢3/4,81 = (U1 — p2)modr and 6 = (u; — p3)mod(2xw/3). If vy =

vzmod 7z then the cosine in (4.14) is equal to —1 and x; = |c; — 3/4 c3|. Hence
the square root in (4.14) is always real. If vi = vsmod2x, then k; = ¢; + 3/4c3,
which is in fact clear from (4.15). O

In this context note that a series expansion for the normalizing constant Gé3)
is provided by (24) in [4]. The precise relationships between the GVMFj and the
GvMy, distributions are straightforward to obtain for the cases iy = 1, k = 1 and
i1 = 1,i3 = 2, k = 2. The assertion that there is an analogue relationship between
GVMF}, and GvMy, densities for 1 < iy < k and k > 3 follows from the fact that
cosk can always be expanded into a sum of multiple angle cosines with multiples
lower than or equal to k. For example, cos® o = (cos 5 4 5 cos 3o + 10 cos o) /16.
Hence, it must always be possible to re-express a GVMF; density (4.1) with 1 <
ir < k in terms of the GvMy density (4.12). However, general formulae for the
GvMy, parameters in terms if the GvMF; parameters seem difficult to establish for
k > 3. So GvMF and GvM distributions are closely related and in fact, Theorem 4.1
and Corollaries 4.1 and 4.2 are the analogues of Theorem 2.1 and Corollaries 2.1
and 2.2 by [4], respectively, of the GvM distribution.

The next results provide characterizations of GvMF distributions in terms of well
known multivariate distributions, Result 4.2 is a characterization in terms of the
multivariate normal distribution. Let us remind that a conditional offset distribution
is the conditional distribution of a random vector given that it has norm equal to one.

Result 4.2. Assume X ~ N (E’ X) in R?, with X positive definite. If I3 # 0, then
X has a GYMF,; conditional offset distribution on S?~!. More precisely,

X|1X] =1 ’\’GVMFPJ,_l(ml,...,mp+1;C1,...,Cp+1;1,2,...,2),

where, given V = (v,,,... ) nonsingular such that ¥~1 = V V7™, we have

%
Vop
my o= [Tl e = 1ZT o myy = el e ¢ =
—||y°j||2/2,f0rj =1,...,p.If p =0, then

XX =1 ~GvMFp(my, ... ,m, 43¢, ..., Cpt152,...,2).

Proof. The conditional random vector X | ||X|| = 1 has density at x € SP~!
proportional to

1
exp ETZ‘_lg—EfZ‘_lg . (4.16)
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Clearly, EZ‘_IK =cimyx. Ifp # 0, then we can define m; and c; as in Result 4.2.
If p = 0, then the term ¢, mx vanishes from the GYMF, 1 density. The three

following statements are equivalent: X is positive definite, X! is positive definite
and, third, there exists V nonsingular such that X~! = VV7. As v, ; denotes the

jthcolumnof V,j =1,..., p,wehave VV" = Zj_l Y Oj With this,

j=1 j=1
p
= ZCJ-H@;'-HE) )
j=1
which terminates the proof. Note that because V' is nonsingular, v,y ....v,, are
linearly independent and the same holds for m,,....m, ;. |

Result 4.3 below extends the characterization of the GvMF distribution given by
Result 4.2 to the situation where the covariance matrix is singular.

Result 4.3. Let X' be a p X p semi-positive definite matrix of rank 7 < p and denote
by A1,...,Ar > 0its non-zero eigenvalues. Let X € R? have the singular normal
density at x € R? given by

r 1
R S (e IR CEy 1) B CA )
where X'~ is a generalized inverse of ¥, i.e., a matrix satisfying ¥ ¥~ Y = X.
Then, for I3 # 0, X has the GYMF,((my,...,m, ic1,...,¢r4151,2,...,2)

conditional offset distribution on S?~!, where the parameters m ;, c;, for j =
1,...,r + 1, have the same form as under Result 4.2, with ¥~ replacing ¥ ~!.
If& = 0, then X has a GYMF, (m,,...,m ;c2,...,¢r+1;2,...,2) conditional
offset distribution on SP~!.

Proof. For A1 = diag(Ay,...,A,) and 7 = (Zol,...,zor), where Yo is the
normed eigenvector of X associatedto A;, j = 1,...,r, wehave X~ = [MAT I}
(see, g, [11, p. 477]). Define V = (vyy.... O,) = NA]Y? Then 3~ =
VVT = Z As the conditional offset density at x € S p 1 is proportional
to

=10 Yo,
Ty — 1T—
expip ¥ z—izﬂz ,

it follows that £" X~ x = ¢ym}x and
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Note that the density (4.17) of X is defined on the translated sub-vector space
{x e R?|N"(x — u) = 0}, where N is a p x (p — r) matrix satisfying N'¥ = 0
and N'N = I,_,. Given ¥ = 'AI'", with A = diag(A1,...,4,,0,...,0) and
I' = (I, I;) with orthogonal and normed columns, X can be expressed as X =
Y + u, where Y ~ N(0, Aq). Hence N can be taken equal to I'>. For further
details refer e.g. to [11, p. 42].

Gatto and Jammalamadaka [5, Characterization 2, p. 348], shows that for the case
p = 2, the conditional offset distribution of a bivariate normal vector is a GvM;
distribution. As noted by Result 4.1 and by the paragraph following it, any GvM;
distribution can be re-expressed as a GvMF, distribution, and hence Result 4.2
generalizes this characterization of the GvM, distribution to dimensions p > 2.
Result 4.2 with u = 0 yields the GvMF,, conditional offset distribution having p
quadratic summands only. In agreement with this, when p = 2, we can see from [5,
Characterization 2], that & = 0 leads to the bimodal von Mises conditional offset
distribution, i.e., to the GVM, density given in (4.12) with k1 = 0.

We can also deduce directly from (4.16) thatif ¥ = 02],foro > 0, and n#0,
then X | || X]|| = 1 ~ vMF(|| ||~ ; 072||||), which is well known.

The next result shows that the GYMF distribution arises also as conditional offset
distribution from the exponential spherical class suggested by [3] as a general-
ization of the multivariate normal distribution. This class consists of multivariate
distributions with densities at x € R? proportional to

exp |~ 1~ W - bt @.18)

where A € R?, W is a p x p positive definite matrix and r > 1.

Result 4.4. Assume X € R? has the exponential spherical density proportional to
4.18) with W = wl, forw > 0,andk =r/2 € {1,2,...}.If A # 0, then X has a
GvMF;, conditional offset distribution on S?~1. More precisely,

X X]| =1~ GvMFi(m,...,m;c1,...,ck;1,...,k),

where m = ||A||~'A and

k

e =2 (M) e @.19)
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forr = 1,...,k. If A = 0, then the conditional offset distribution of X is the
isotropic one.

Proof. The conditional random vector X | ||X]|| = 1 has density at x € SP~!
proportional to

wk
exp § =[x = 1) (x - )
The above exponent can be expanded as

r k

w k T _N\F Ty \k—r
—_ -2 1+
2k,=o(r)( Ax) (1 + A7,

which is, up to an additive constant and for A # 0, equal to Zlle cr(m™x)", with
m and ¢, as given above, forr = 1,... k. O

From Result 4.1, the GvMF; distribution can be reparametrized in the GvM3
form when p = 2 and thus Result 4.4 extends in some way the characterization
of the GvM, distribution in terms of the bivariate normal distribution given in [5,
Characterization 2]. The case k = 3 is given precisely in Result 4.5 below.

Result 4.5. Assume X € R? has a density proportional to (4.18) with W = w1, for
w > 0and k = r/2 = 3. Then

arg{ X} [ ||X]|| = 1 ~ GVvM3 (1, ph2, U3 K1, k2,K3),

where for c;, ¢p and c3 given by (4.19), u; is given by (4.13) and k; by (4.14) with
vy = v3 = arg{A}, no = u3 = arg{r}, ko = ¢3/2 and k3 = c3/4.
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Chapter 5
A New Nonparametric Test of Symmetry

Kaushik Ghosh

Abstract We present a new nonparametric test for symmetry about an unknown
location and investigate its large sample properties. Asymptotic normality of the test
statistic is established and an estimator of the asymptotic variance is also presented.
Results of a simulation study and data analysis are also presented.

5.1 Introduction

In many practical situations, it is important to determine whether the underlying
population has a symmetric distribution. For example, if one is interested in esti-
mating the measure of location, having a skewed distribution would give rise to
consideration of more than one such measure. In the case of a paired sample prob-
lem with “treatment” and “control”, the hypothesis of “no treatment effect” implies
that the paired difference Z = X — Y is symmetric about 0. Many robust statisti-
cal methods (see [35]) depend on the assumption of symmetry. In case symmetry
is not valid, one would need to determine a symmetrizing transformation before
applying the statistical procedures. Koziol [40] mentions using a test for symme-
try as a screening procedure before applying the modulus family of transformations
introduced by [36] for bringing data to be closer to normality.

The problem of testing univariate symmetry has received attention in the lit-
erature for quite some time. Past work in this area can be broadly classified
into two groups — one where the center of symmetry is known (without loss
of generality, this is then taken to be 0) and the other case is where the cen-
ter of symmetry is completely unknown. A common class of nonparametric tests
in the first category is the weighted sign test, which has been studied in detail
by many authors, such as [29, 30, 37, 59]. Special cases of the former include

K. Ghosh
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the popular sign test and the signed-rank test. Other examples include works by
[14,20,47,53,56]. Aki [3] proposed a new test of symmetry which generalized the
results of [14] and [53]. Ahmad [1] proposed asymptotically normal estimates of
symmetry statistic by [53]. Reynolds [51] and Rosenstein [52] developed a sequen-
tial signed-rank test for symmetry about a known value. Further work have been
done by [6,8,9,17,21,31-33,39,41,42]. Some of the recent work in this area have
been by [2, 15,45, 46, 58].

The case of unknown center of symmetry is a more realistic and practical prob-
lem. One of the earliest articles in this area is by [34] where the authors use the
difference of the mean and median as a measure of skewness. One group of tests is
obtained by applying the tests with known center, where the center is estimated from
the data. Gastwirth [26] proposed the modified sign test, whereby the unknown point
of symmetry is estimated by the sample mean and a sign test is developed for sym-
metry around this point. Other articles in this category include those by [10,21,28].
Boos [13] propose a test for symmetry based on the Hodges—Lehman estimator of
location. Davis and Quade [19] and Randles [50] propose asymptotically distribu-
tion free tests of symmetry. Antille and Kersting [5], Antille et al. [6] and Finch [25]
propose tests based on symmetric differences of spacings. Csorgd and Heathcote
[18] and Koutrouvelis [38] use the empirical characteristic function to develop tests
for symmetry about an unknown location, that do not require estimation of the
unknown location. Koziol [40] propose using rank-based tests of symmetry with
known center where the center is estimated using asymptotically efficient methods.
Eubank et al. [23] also propose a class of rank-based tests of symmetry. Schuster and
Barker [54] proposed a symmetric bootstrap procedure for testing symmetry about
unknown center, which was later examined in further detail by [7] and extended to
the dependent case by [48]. Cabilio and Masaro [16] propose a test of symmetry
about an unknown location based on deviation of the sample mean from the sam-
ple median, similar to the lines of [34]. Some of the recent work in this area are
[22,43,44].

In this article, we develop a new test of symmetry for univariate distributions
when the center of symmetry is unknown. Our test is based on a new characteriza-
tion of symmetry. This article proceeds as follows. In Sect. 5.2, we propose the new
estimator and investigate its properties. In Sect. 5.3, we provide results of simulation
studies in support of the theoretical properties. In Sect. 5.4, we provide a real-life
example of use of the proposed statistic. Finally, we end with concluding remarks
in Sect. 5.5. The proofs of results presented here are presented in the Appendix.

5.2 Theory

Suppose X7y, ..., X, is a set of observations (not necessarily i.i.d.) from an under-
lying continuous distribution with cdf F' and corresponding pdf f. We want to test
for symmetry of f about an unknown point u (say). For any p € (0, 1), consider
the function
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F ' (p)+F'(1 —p))_

g(p)=F( 7

Note that g(p) < %iff is left skewed, g(p) > %iff isright skewed and g(p) = %

is f is symmetric. Also note that g(p) = g(1 — p). Consequently, we propose the
following quantity to characterize the symmetry of f:

1/2
r=t(F) = /0 ¢(p)dp. 5.1)

It follows from prior remarks that t = i for symmetric, t > i for right skewed and
T < % for left skewed distributions.
Suppose Fisan empirical-process based estimate of F', such that

N 2 (5.2)

where WF is a Gaussian process with covariance kernel Kfr(-,-). We propose
estimating t using the quantity

1/2 —1 r—101 _
%:/ F(F P+ F 0 p))dp. (5.3)
0

2

Our goal is now to estimate the asymptotic distribution of 7. Consider the map
¢ : D[0, 1] — DJ0, 1] given by

(5.4)

F'(p)+F (1 —p))
5 .

¢(F)(p) = F (
We first present an important result.

Theorem 5.1. Suppose F is a distribution with pdf f and F be an empirical-
process based estimate of F satisfying (5.2), where WF is a Gaussian process with
covariance kernel K (-, -). Suppose we define the functional G = ¢(F) and G be
its empirical counterpart, where ¢ is given by (5.4). We then have

V(G -GS wg.

where Wg is a Gaussian process with covariance kernel Kg(-,) given by

Ko(s.1) = Kr (F_‘(S) +§—1(1 -5 F0) +§_1(1 _t))

1 (F Y s)+F'(1-ys) 1 _ F ')+ F~1(1-1)
-/ 2 ) 7 e (o 2 )
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ﬁ ( S, _1“”;““(1—0)}

_§f( 1<t)+2F 1(1—t)){fF11(t) ( o _I(S)+2F—1(1_s))
+ﬁm (F—1(1_,), 1(s)+§—1(1_s))}
+%f(F—‘(s)+§“(1—s))f(F—‘(t)+§—1(1_t))

X{ K (F@. F()

FF1(s) FF1(1)
! F(F1(1—5), F71(1))

e

1 - -1
+fF—1(s)fF—1(1—t)KF (F~'(s). F~'(1—=1))
1

T =Y =T

F(F'(1—s), F7'(1— t))} .

Corollary 5.1. Suppose X1...., Xy, areiid. F, which is symmetric about u and
F is the corresponding empirical distribution function. We then have

t }+1 )
JF~ 1() fE1@) 2fF () fF71()

Theorem 5.2. Suppose X1, ..., X, are observations from a distribution F. Let T
and T be as defined earlier. We then have

Ka (s, t)—— f( ) (s AT).

Vi —1) 5 N, 0?)

where

1/2 p1/2
w? = / K (s, t)dsdt.
0 0

Corollary 5.2. Suppose X1,..., X, are i.i.d. F which is symmetric about ji. Fur-
ther, assume that F has finite second moment. We then have

Jn (% - %) LN, 0?),

where |
o = E——f(M)E(IX b + f (WEX — ). (5.5)
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Table 5.1 Asymptotic variance of the symmetry statistic for some special symmetric distributions

Distribution w?
Uniform ‘% = 0.0208
Normal ”1—2 = 0.0227
Laplace + =0.0625
2
A ret s
t(v>2) i { r(g) } o= De=D)
2
1 m—+1 r(2m)
Beta(m’ m) 16 16"m(2m—-1) {Fz(m)}
— 2
Logistic % = 0.0273

Note that the expression for w? is invariant with respect to location and scale

changes. Also note the similarity of the asymptotic variance formula to that of the
asymptotic variance of Sk defined in [16].

Table 5.1 gives the asymptotic variance of the symmetry statistic for some special
distributions. It is interesting to note that the asymptotic variance for the case of
samples from ¢, distribution decreases with v and — == —2 a5 v — oo. Similarly,
for samples from the Beta(m, m) distribution, the asymptotic variance — ’lrﬁ—n as

m — oo and — E as m — 0. The minimum is attained at m = 1.66529.

5.2.1 Estimation of the Asymptotic Variance

The asymptotic variance w? of the 7 statistic given in (5.5) depends on unknown
population quantities, which need to be consistently estimated. From [57] and [12],
we see that the variance of 1 37 | |X; — X| is of bounded order O(n™") and
ELST [ 1Xi — X| = E|X — pl{l + O(n™/2)}. Hence, 1 3°7_ |X; — X is
a consistent estimator of E|X — u|. Consequently, we can consistently estimate w
along the lines of [16] using

o* = ———f() ZIX Al+ 7 f(M)Sz (5.6)

1—1

where S? = ﬁ (X — X)? is the sample variance. Here [ is the sample
median and f (f0) is the kernel density estimator of f (i) obtained using

1 & x—X;
nhnEK( hy, )

where K(-) is the Epanechnikov kernel

fx)=

3 1
K(t) = { 7z (1= 517 for || < V/5
0

otherwise,
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and the bandwidth #,, is chosen to be
h, = 1.06 min(std. dev, IQR/1.34)n"'/3

(see [55, pp. 43—47] for more on these choices).
Note that for a given random sample X1, X», ..., X, we can write

%[#<X5m> +#(X5%) +...+#(X < M@)] if n is even

Ll (x < 2oti) gy (x < dotfemn) 4y g (X<Xon )] ifnisodd

Hence, a 2-sided test for symmetry at level of significance o can be given as:

reject Hy if Z = \/n

s 1
Iz
w

> Za/2-

5.3 Simulation Studies

We conducted a simulation study based on samples generated from a generalized
lambda distribution (GLD) considered in [42, 58]. The quantile function of the
generalized lambda distribution is given by (see [49])

uts — 1- u)M

x=Fl'u) =1+ P ,

0<uc<l.

Here A7 is the location parameter of the distribution, A, the scale parameter and
A3, A4 determine the skewness, with symmetry when A3 = A4.

Data were generated from 14 members of the GLD family. Foreach (A1, A2, A3, A4)
combination, we considered 4 sample sizes (n = 30, 50, 100 and 500). For each
n, we generated a random sample of size n and performed four different tests of
symmetry about an unknown location: (i) the proposed test (denoted by 7), (ii) the
test proposed by [43] (denoted by T'), (iii) the test proposed by [44] (denoted by y1)
and (iv) the test proposed by [16] (denoted by C).

Note that each of the 4 test statistics are asymptotically normal whose asymp-
totic variances depend on the underlying distribution. In practice, one may decide to
estimate this asymptotic variance along the lines of Sect.5.2.1 to make these tests
completely distribution free. However, a brief simulation study (not presented here)
indicated that the resulting tests have poor behavior, especially for small samples.
This is possibly due to the slow convergence of the estimator of the asymptotic
variance and/or due to slow convergence to normality.

Consequently, we decided to follow the methodology used by [16, 43, 44],
whereby the critical values of the 4 tests are first empirically estimated based on 10°
simulations from a normal distribution. The resulting critical values are then used to
study the power of the tests, with each power calculation based on 10° simulations.
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Table 5.2 Estimated type I error probabilities of the various tests of symmetry when data are
simulated from some symmetric members of the GLD family. Critical values are based on normal

reference distribution. Results are based on 10° simulations, with nominal & = 0.05

Test P(type I error)
n =30 n =50 n = 100 n = 500
Case 1: A = (0,2,1,1)
T 0.0443 0.0495 0.0437 0.0411
T 0.0940 0.1029 0.1006 0.1064
Vi 0.1151 0.1271 0.1308 0.1376
C 0.1173 0.1295 0.1314 0.1376
Case 2: 1 = (0,0.19745,0.134915, 0.134915)
T 0.0539 0.0498 0.0507 0.0476
T 0.0531 0.0523 0.0518 0.0501
Y1 0.0524 0.0491 0.0515 0.0496
C 0.0541 0.0505 0.0521 0.0496
Case 3: A = (0, —1,—0.08, —0.08)
T 0.0928 0.0930 0.0938 0.0942
T 0.0609 0.0625 0.0635 0.0588
Y1 0.0409 0.0446 0.0433 0.0382
C 0.0421 0.0457 0.0439 0.0381
Case 4: A = (0,—0.397912, —0.16, —0.16)
T 0.1106 0.1229 0.1178 0.1344
T 0.0806 0.0827 0.0769 0.0773
Vi 0.0459 0.0446 0.0444 0.0439
C 0.0476 0.0463 0.0449 0.0438
Case 5: A = (0,—1,—0.24, —0.24)
T 0.1416 1475 0.1585 0.1820
T 0.1016 1069 0.1061 0.1074
Y1 0.0457 0.0475 0.0454 0.0491
C 0.0478 0.0482 0.0461 0.0491
Case 6: A = (0,—5.11256, —1, —1)
T 0.4300 0.4771 0.5413 0.6621
T 0.5364 0.5908 0.6663 0.7983
Y1 0.0945 0.0864 0.0907 0.0991
C 0.0987 0.0895 0.0912 0.0989
Table 5.2 gives the results for the symmetric cases (A; = A,). We see from

Table 5.2 that for case 1 (uniform), the only recommended test is 7, since it is the
only one maintaining the type I error level. For cases 3, 4 and 5, tests 7 or T should

not be used — instead one should preferably use y;. For case 6, none of the four
tests are recommended. Finally, note that all the tests more or less maintain their

significance levels for case 2. This is expected, since case 2 gives rise to a normal-

like distribution.

Table 5.3 gives the results for the asymmetric cases. The density curves of the dis-
tributions are sketched in [42] and denoted there as case 2 to case 9. From Table 5.3,
we see that for case 11, all the tests have very low power, even for quite large sample
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Table 5.3 Estimated power of the various tests of symmetry when data are simulated from some
asymmetric members of the GLD family. Critical values are based on normal reference distribution.

Results are based on 10° simulations, with nominal « = 0.05

Test Power

n =30 n =150 n = 100 n = 500
Case 7: A = (0,1,1.4,0.25)
T 0.1872 0.2759 0.4627 0.9884
T 0.2692 0.3756 0.5668 0.9908
Y1 0.2808 0.4002 0.5993 0.9933
C 0.2851 0.4041 0.5994 0.9933
Case 8: A = (0,1,0.00007,0.1)
T 0.6048 0.8141 0.9812 1.0000
T 0.6563 0.8388 0.9815 1.0000
Y1 0.5972 0.8111 0.9787 1.0000
C 0.6032 0.8145 0.9788 1.0000
Case 9: A = (3.586508, 0.04306, 0.025213, 0.094029)
T 0.2482 0.3850 0.6368 0.9993
T 0.2375 0.3533 0.5952 0.9985
Y1 0.2019 0.3201 0.5690 0.9984
C 0.2071 0.3256 0.5696 0.9984
Case 10: A = (0,—1,—0.0075, —0.03)
T 0.3981 0.5979 0.8678 1.0000
T 0.3793 0.5748 0.8424 1.0000
Y1 0.3035 0.5001 0.8058 1.0000
C 0.3107 0.5063 0.8064 1.0000
Case 11: = (—0.116734,0.351663, —0.13, —0.16)
T 0.1350 0.1489 0.2021 0.5145
T 0.0995 0.1076 0.1339 0.4132
Y1 0.0565 0.0636 0.0879 0.3290
C 0.0587 0.0648 0.0885 0.3285
Case 12: A = (0,—1,—0.1,—0.18)
T 0.2628 0.3813 0.6058 0.9974
T 0.2224 0.3279 0.5345 0.9927
Y1 0.1309 0.2217 0.4249 0.9874
C 0.1360 0.2259 0.4255 0.9873
Case 13: A = (0,= —1,—0.001, —0.13)
T 0.7935 0.9490 0.9989 1.0000
T 0.8398 0.9624 0.9986 1.0000
Y1 0.7545 0.9370 0.9981 1.0000
C 0.7608 0.9390 0.9981 1.0000
Case 14: . = (0,—1,—0.0001, —0.17)
T 0.8260 0.9616 0.9998 1.0000
T 0.8724 0.9715 0.9999 1.0000
Y1 0.7908 0.9491 0.9997 1.0000
C 0.7960 0.9502 0.9997 1.0000
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sizes. For cases 9, 10, 11 and 12, the test T seems to be slightly preferable over the
other choices. Note that in all these cases, the distribution is close to symmetric and
is thus hard to distinguish. In none of the 8 cases, the test y; seems to be preferable.

5.4 An Example

We use the data from [20] to illustrate our proposed method. Table II of [20] (repro-
duced in Table 5.4) gives survival times (in days) of 72 guinea pigs that received a
dose of tubercle bacilli (see [11] for more details on the data). A plot of the kernel
density estimate of the data is given in Fig.5.1.

Table 5.4 Survival times (in days) of guinea pigs. Data reproduced from [20]

10 33 44 56 59 72 74 77 92 93
96 100 100 102 105 107 107 108 108 108
109 112 113 115 116 120 121 122 122 124
130 134 136 139 144 146 153 159 160 163
163 168 171 172 176 183 195 196 197 202
213 215 216 222 230 231 240 245 251 253
254 254 278 293 327 342 347 351 402 432
458 555

Estimated density of survival times of 72 guinea pigs

Density
0.003 0.004 0.005
1 1 1

0.002
1

0.001
1

0.000
1
|
\

T T T
0 200 400 600
Days

Fig. 5.1 Kernel density estimate of the guinea pig survival times
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Application of the proposed test resulted in a test statistic value of Z =
1.597538, which corresponds to a p-value of 0.0550 against the alternative hypoth-
esis of positive skewness. Thus, the data is close to significantly positively skewed
at 5% level.

5.5 Conclusion

We have proposed a new test of univariate symmetry about an unknown loca-
tion. From simulation studies, the test is seen to possess improved power over
other choices when the underlying distribution is close to symmetric. The results
presented here are based on estimating the critical values assuming a normal distri-
bution. Preliminary simulations have shown that the asymptotically distribution-free
version (obtained by estimating the asymptotic variance) has poor performance,
especially for small sample sizes. We believe that this is due to the slow conver-
gence to the limiting distribution. Methods to speed up convergence need further
investigation.

Although we have presented the expression for the asymptotic variance only for
the case of i.i.d sample, the same principle can be used for cases where one has
ranked set samples or its generalizations (see [27]). The case of testing symme-
try based on such generalized sampling schemes is currently ongoing and will be
presented in a later article.

Appendix

Lemma 5.1. Consider the map ¢ : D[0, 1] — DI0, 1] given by

sy = F (L2 AED),

2

Let ¢ (h) be the Hadamard derivative of ¢ (F) in the direction of h. Then, we have

-1 1 _ 1 1 B
e e 2 R

y [h(F_‘(p)) n h(F~'(1 —p))}
SFEp)  fFETA=p)]

Proof of Lemma 5.1: The Hadamard derivative of ¢(F) in the direction of 4 is
given by
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¢ (F +th)(p) — (F)(p)
t

b (1) (p) = lim

= lim ~[$(F + 10)(p) ~ $(F)(p)]

_ }E‘},% [(F i) ((F + th)~(p) +§F + )71 — p))
_F(F“(p)+f“(1—p))]

1 (F 4+ th)y Y (p) + (F +th)"'(1 — p)
Z}%?[F( 2 )
e ((f + th) "' (p) +§F + th)~' (1 — p))

_F(F“<p)+f—1(1—p>)'

, 1[ ((F+th)_1(p)+(l_7+th)_1(1—p))
=lim-|F

t—0 2

_F(F_I(P)"’;_l(l—P))_

+h((F+th)_1(p)—i—;F—}-th)_l(l—p)).

Letu = F- Y (phu+v = (F+th Y (pha=F'1l-p.a+b=(F+
thy™'(1 — p). Then, Fu) = p = Fu+v) +th(u+v)and Fla) = 1—p =
F(a+ b) +th(a + b). Note thatas t — 0, u — 0 and v — 0. Hence,

N ((F +th)H(p) + (F +h)~'(1 —p)) _ (u+v+a +b)
2 2

u-+a
—>h( 5 )
_ h(F‘l(p)JrF‘l(l—p))
5 )

(5.7)

Also,

1[F((F +th~'(p) + (F +th)7'(1 —p)) _F(F_l(p) +F71 —P))}
t 2 2

u+ v+b u+
F(Ta+T)_F(Ta) b
E z

=T} x T5 say.
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Ast — 0, we have

o f (u _; a)
-1 —101 _
_ f(F (p)+;F a p)). (5.8)
Also,ast — 0,
p Y +b
Y
1 vh(u +v) bh(a + b)
T2 |:F(u) —F(u+v)  F(a)— F(a+ b)}
N l (—h(u) n —h(a))
2\ fw)  fla)
_ —L[hF(p) | h(F7'Q —P))]
-2 [f(F“(p)) N GErE G2
Combining (5.7), (5.8) and (5.9), we get the result. O

Proof of Theorem 5.1: From (5.2) and the functional delta method (see [4,24]), we
have

Ji@(F) = $(F)) S ¢ (Wr) (5.10)
where
-1 —101 _
$r (W) = wr (L)
Ly (F‘l(p) +F7'1 —p))
2 2
§ [WF(F—‘(p» LW —p))}
fF=Y(p)) fF1d=p) |
The result follows from the above. O
Proof of Theorem 5.2:
1/2 . 1/2 d 1/2 p1/2
ﬁ( ¢(F)(p)dp —/ ¢(F)(p)dp) —>/ Kg (s, t)dsdt.
0 0 0 0

Proof of Corollary 5.2: Note that
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1/2 p1/2 s 1 [#
/0 /0 fF_—l(s)dsdt = 5/_oo(u—t)f(t)dt-

Also,
1/2 p1/2 SAL m
dsdt = / (w =12 f(@t)dr.
/o o JFU)SF() —o0
Combining, we get the result. O
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Chapter 6

A Semiparametric Bayesian Method
of Clustering Genes Using Time-Series
of Expression Profiles

Arvind K. Jammalamadaka and Kaushik Ghosh

Abstract An increasing number of microarray experiments look at expression lev-
els of genes over the course of several points in time. In this article, we present two
models for clustering such time series of expression profiles. We use nonparametric
Bayesian methods which make the models robust to misspecifications and provide
a natural framework for clustering of the genes through the use of Dirichlet pro-
cess priors. Unlike other clustering techniques, the resulting number of clusters is
completely data driven. We demonstrate the effectiveness of our methodology using
simulation studies with artificial data as well as through an application to a real
data set.

6.1 Introduction

Rapid advances in genome-sequencing technology has made it possible to collect
data simultaneously on expression-levels of all genes of an organism relatively eas-
ily than it has been ever before. Usually, the number of genes is in the thousands
and the data might be very high-dimensional, thus requiring specialized statistical
tools for analysis. One of the natural questions that scientists have been pondering
over is how to use such expression data to cluster genes into functionally related
groups. The aim is thus to identify genes with similar expression patterns, which
can further lead to improved understanding of cell functioning — possibly opening
way for better treatment of diseases.

The expression data can be in the form of a single snapshot at a point of time or
can be in the form of multiple measurements of the genes, taken at several points
in time. The latter gives rise to time series of expression profiles and is naturally
more informative in clustering genes than if one were to use single time snapshots.
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Time series experiments are becoming increasingly common (see [2,8-10,12,13,15]
for some recent examples of work in this area) with the improvements in high-
throughput data collection techniques such as microarrays.

Analyzing such expression data present several special challenges, since one
needs to account for the correlation among several genes at a timepoint as well as
correlation among measurements on the same gene at different timepoints. Several
of the past methods dealing with such data have assumed that the expression profile
for each gene is a multivariate vector where the profile vector, given the parameters,
is assumed to be multivariate normal with independent components (see, e.g., [8—
10]). While such simpler models provide a first-cut, they fail to take into account the
dependence of the measurements on a gene at various timepoints and are thus not
realistic. In this paper, we in contrast, allow for potentially rich correlation struc-
tures between timepoints. Such dependence between expression values for the same
gene, is quite natural in our opinion, and is the main contribution of this paper.

This paper is organized as follows. In Sect. 6.2, we present two proposed models
for the time series of expression data. In Sect. 6.3, we discuss the techniques used to
estimate the model parameters. To demonstrate the ability of the proposed models
to correctly identify the underlying clusters, we present results of a short simulation
study in Sect. 6.4. In Sect. 6.5, we illustrate this method by an application to the
well-studied yeast cell-cycle data [14]. Finally, we present concluding remarks in
Sect. 6.6.

6.2 Model Description

Suppose that a microarray experiment consists of N genes. Each gene is repeat-
edly measured over time on p different occasions, resulting in a p-dimensional
measurement vector y; = (y,-l,...,y,-p)’ forgenei (i = 1,...,N). Let u; be
the mean vector of y; and X; be the corresponding covariance matrix. In what
follows, we present two different models for the joint distribution of the measure-
ment vectors y;. The models differ in how the variance-covariance matrix X'; is
represented.

6.2.1 Heteroscedastic Model

In this model, we assume that the variance-covariance matrix for the measurements
on gene i is givenby X; = UI-ZH (pi). Here 01-2 and p; are gene-specific variance and
one-step correlation parameters respectively and H(-) is a matrix-variate function
that is common to all genes. The matrix H(-) provides the dependence structure
among the measurements at various timepoints on a particular gene and is assumed
to be completely known.
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Two possible forms of the H(-) matrix are

H(p) = (0" 1)) pxp

and
H(p) = ((Li=j3 + pLii#53) pxp-

The first form assumes that the correlation between measurements at any two
timepoints decays exponentially as a function of the distance between the two time-
points, so points further away are less correlated with one another. The second form
assumes that the correlation remains the same between any two points of time.

We assume that conditional on the mean vector, the common variance and
one-step correlation, gene expression profiles for various genes are independent
multivariate normal random vectors. That is,

indep

yil(rioofs pi) "~ Np(uis o7 H(pi)),  i=1,....N.

Let 0; = (u;, 07, p;) be the vector of parameters used to describe the joint
distribution of the expression profile of ith gene. We will say two genes i and j
cluster together if their corresponding parameter vectors #; and 6 ; are identical. To
this end, we assume that 6; |G % G and G|(M,Gy) ~ D(M, Gy). Here D(M, Gy)
denotes a Dirichlet process [5] with base measure G and concentration M .

The reasons for such a choice of prior distribution are 2-fold. First, using a
Dirichlet process makes the distribution of the parameter vectors flexible and robust
to mis-specifications. Second, due to the natural clustering property of samples
from a distribution with a Dirichlet process prior [1], there is inherent data-driven
clustering among the model parameters.

We assume that under the baseline prior Go, (1;, 67, p;) are independent with
multivariate normal, inverted-gamma and beta distributions, respectively. In other
words,

Go = Np(po, Xo) x IG(ag., bs) x Beta(a,, by)

where

Yo = diag(og,, . . . ,ng)
with

02,02, X 1G(ao, bo)
and

o = (o1, .- wMOp)T-
We further assume

M ~ Gamma(ap,bpy).

All the hyperparameters aq,bg,a0,bo,ap,bp, am.bm, o1, ..., lop are
assumed known and in practice, are chosen to make the priors as noninformative
as possible. Due to lack of closed form expressions for the posterior distribution
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and easy availability of univariate conditionals, we will use a Markov chain Monte
Carlo approach to estimate the model parameters. The natural clustering property of
the Dirichlet Process will lead to clustering of the values of ;.

6.2.2 Homoscedastic Model

In this case, we assume the variance of measurements is same across all genes and
across all timepoints. Hence, the clustering of genes is achieved based on the gene-
specific mean vector and one-step correlation. Thus, we have

inde, .
yil(u;. 02 pi) "~" Np(u;.02H(p;)). i=1,....N,

where N = number of genes,

H(p) = (0" pxp,

or
H(p) = ((Igi=jy + pli#j3) pxp-
is the correlation matrix, and p = number of observations per gene.
We further assume:

iid
(ki,pi) =06 ~ G,

G ~ DP(M, Gy),

Go = Np(pg, Xo) x Beta(ap, bp).

Yo =diag(o}, ,olz,)pxp,
0r.....0, “1G(a*, b*),

Ro = (o1, - -, Hop)'s

M ~ Gamma(ap,bar),

0% ~ IG(ag, bs).

Note that this model differs from the heteroscedastic model by the fact that all
genes are assumed to have the same variability in their pointwise measurements and
cannot be obtained as a special case of the former model.

6.3 Model Fitting and Estimation

Due to lack of closed-form expressions of the posterior distributions, model fitting
is done using Markov chain Monte Carlo procedures. In particular, since univariate
conditionals of the model parameters are easily obtained, we use the Gibbs sampler.
Details of the implementation of the Gibbs sampler are outlined below.
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6.3.1 Heteroscedastic Model

We can update the model parameters according the following scheme:

-1

N 1 1 N
2 2
00j|"""1G ao+7,|:a+§;1(ﬂij_ﬂoj):| s

j=1...,p.

2. Due to the clustering property of samples generated from Dirichlet process,
not all values of 6; are distinct. In any iteration, let there be k distinct values,
0F,....0;. Let¢ = (c1,...,cn) be the configuration vector where ¢; = j if
and only if 6; = 9}‘, Jj = 1,...,k. Let n; be the number of occurrences of 9}‘.
The configuration vector is updated every iteration using algorithms of [11].

3. Update the distinct values 9;-‘ as follows (j = 1,...,k)

2 2
fasor o5l o [T fOilisof .05 f(uhlio. Zo)

ici=j
2
x f(0} lac.bs) f(pjlap. by).
Consequently, it is easy to show that

@ |-+ ~ NGrj, I}f) where

-1

*2 -1
re=| (2 mpy) +5
i = " Pj 0

J
and
o - 1
=T} (#H(p}f)) — 2 v+ e
(b) J T ;=
-1
2 pn; 1 1 _
o |-~ 1G aa+T’,[b—+5 > i —uHTH(Y) I(Yi_ll-j):| .
o . .
ici=j
(c)
1 —L‘*zZ,-;Ci_,(yi—ujf)TH(pj)“(yi—ujf)}
fojl) X———¢ 7
|H(Pj)| 2

x (P (1= pp)tet. 6.1)
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The components of 9;‘ can be updated one at a time using the above results.
Updating of ,o}‘ using (6.1) can be done using Adaptive Rejection Sampling
(ARS) or Adaptive Rejection Metropolis Sampling (ARMS) (see [6, 7]).

4. M is updated using a mixture of Gammas as outlined in [4].

6.3.2 Homoscedastic Model

The parameter updates are done according to the following scheme:

1. Update the pointwise variance using

np | 1 1Y -
2 s N1y, — s

o]~ IG a0+7’|:z+§§(y' wi)H(pi)~" (yi Hz)i|

2. Update the entries of the baseline covariance matrix Xy using

-1

N |1 1g
2 2
oyl ~1G a*+?,[ﬁ+§;1(uu—uw)} :

j=1...,p.

3. As in the heteroscedastic model, let there be k distinct values, 67, ..., 9,:‘ and
let ¢ = (cq,-..,cn) be the configuration vector where ¢; = j if and only if
0, = (9;‘, Jj = 1.... k. Let n; be the number of occurrences of (9;‘. We update
the configuration vector using algorithms of [11].

4. Update the distinct values as follows:

wilo~ Np(u3. I},

o? - !
ry = [(n—j_H(p») ' 2&‘}

where

and

o? 1 _
wi=Tr; (7H(pj)) — > i+ 55 e
J ici=j
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1

f(P;f| ) X ;.
\H(p|

% e—[z(%z Yitej=i Wi —Mj)TH(Pj)_I(J’i—Mj)](pj)ao—l(l _ pj)bp—l_
Use ARS or ARMS to do the last update.
5. Update M using a mixture of Gammas, along the lines of [4].

6.4 A Simulation Study

Performance of both the heteroscedastic and homoscedastic models were evaluated
using a simulation study involving a synthetic data set. This data consists of expres-
sionon N = 60 genes measured at p = 10 equally-spaced timepoints, without loss
of generality which are takentobe t; = 1,...,f19 = 10. The 60 genes are divided
into 3 groups, each consisting of 20 genes. The data were generated according to
the following model
Yij = ¥i(t)) + ey
where
Vi(t) = 8io + 8irt + Bir(t —t1)4 + -+ + Bip(t — 1)+

is the mean trajectory of the ith group and e;; is the random error in measuring the
expression level for gene i at timepoint ;. The parameters for the mean trajectories
for the three groups are given in Table 6.1. The errors for each group are assumed to
be normal with zero mean and variances given by 0.25, 1.0 and 0.04 respectively. In
addition, we also assumed that there is a correlation of p = 0.3 between time-points.

Once we generated the data, the Gibbs sampler was used to sample from the pos-
terior distribution for the fitted models. Both the homoscedastic and heteroscedastic
models were fitted to the simulated data. A burn-in of 10,000 iterations was found to
be enough to reach steady-state. An additional 10,000 iterations were used for the

Table 6.1 Synthetic data trajectory parameters

Group 1 Group 2 Group 3
8io 0 0 —1.0
8i1 0.5 0.5 0.2
Bii 0 0 0
Bir 0 0 0
Bis 0 0 0
Bia 0 0 0
Bis 0 —1.5 0
Bio 0 0 0
Bi 0 0 0
Bis 0 0 0
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Fig. 6.1 Heatmap of clusters of the synthetic data based on the average incidence matrix from
the heteroscedastic model. Higher intensity (in deeper red) denotes more likelihood of clustering
together, while lower intensity (in lighter yellow) denotes less likelihood of clustering. Note that
the three original groups of 20 units each are fully recovered (denoted by the three large squares
on the diagonal)

purposes of posterior inference. For each iteration /, we obtained a configuration
vector ¢ = (cfl), . ,cl(\l,)) of the cluster-structure of the genes. This gave rise to

an incidence matrix Qj(\l,)x N> Where o0 = ((ql.(,l]).)) and ql(l]) = lif cl.(l) = cﬁl), 0

otherwise. The average incidence matrix Q = % ;}1{’ 00 0" was then computed.
The heatmap routine in R was used on 1 — Q to generate a heatmap of the
clustering of the genes based on this average incidence matrix. The result for the
heteroscedastic model is shown in Fig. 6.1.

We note that these models generally seem to perform very similarly on the test
data. While both of them are able to isolate members of the same original group into
clusters, the clustering is not perfect in all the iterations.
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6.5 Application to Spellman Data

The proposed models were next applied to a standard subset of the well-known data
on time series of gene expression of yeast cells due to [14]. Details on the data and
how to access it can be obtained from http://genome-www.stanford.edu/cellcycle.
The subset consists of observations on N = 798 genes measured at p = 18 time-
points. Both the homoscedastic and the heteroscedastic models were fit to the data.
A burn-in of 10,000 iterations stabilized the sampler. After that, an additional 5,000
iterations were used to estimate the model parameters. Each iteration produced a
configuration structure of the genes that cluster together, giving rise to an incidence
matrix. The average incidence matrix was used to ascertain the “average cluster
structure” of the genes using the method of [3] or through heatmaps as described
earlier. The resulting heatmap for the heteroscedastic model is provided in Fig. 6.2.

Gene #

Gene #

Fig. 6.2 Heatmap of clusters of the 798 genes in the Spellman data based on the average incidence
matrix for the heteroscedastic model. Higher intensity of red denotes more likelihood of clustering
together
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Fig. 6.3 Histogram of number of clusters generated using the heteroscedastic model on the
Spellman data
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Fig. 6.4 Mean curves of top 5 clusters found by applying the heteroscedastic model to the
Spellman data
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The median number of clusters using the homoscedastic model is 5 with a 95%
credible interval being (1, 13) and the corresponding figures for the heteroscedastic
model were 7 and (2, 14) respectively. The histogram of the number of clusters in
the heteroscedastic model is given in Fig. 6.3. All the above models were coded in
C, and the code can be obtained from the authors upon request.

A plot of the mean expression profiles for the top 5 clusters corresponding to
the heatmap and the resulting dendrogram for the heteroscedastic model is given in
Fig. 6.4. To get a rough measure of cluster separation, we consider a rather crude
measure of distance achieved by each of the (cluster-specific) average trajectories
from the overall mean trajectory y, as measured by the sum across the 5 clusters and
the 18 timepoints using

5 18
DO i —=)>
i=1t=1
This measure has a value of 39.68 for the heteroscedastic model and 40.13 for the
homoscedastic model. Hence, in terms of the measure of separation, of the clusters
the two models are quite close.

As a check of how robust our choice of prior parameters on M are, we changed
these to yield an expected number of clusters that is closer to 5. The resulting mean
curves for the top 5 clusters are nearly indistinguishable from those obtained previ-
ously. This assures us that the hyperparameters do not play a very significant role,
which is often a point of concern in Bayes methods.

6.6 Conclusion

We have provided a semiparametric Bayesian approach to clustering genes based
on their expression profiles, which we assume have certain correlation structure.
This method is easily implemented using standard computer languages and produces
results in a reasonable period of time (it took about 2 hours to cluster the 798 genes
on an Apple MacBook Pro with 2.6 GHz Intel Core 2 Duo and 4 GB of RAM). One
can easily add additional steps to the Gibbs sampler to make the model more robust
to mis-specifications (e.g., using a multivariate ¢ instead of multivariate normal).
The model can also be used to handle missing data by replacing them with their
imputed values or as an additional MCMC step.
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Chapter 7

On Implementation of the Markov Chain
Monte Carlo Stochastic

Approximation Algorithm

Yihua Jiang, Peter Karcher, and Yuedong Wang

Abstract The Markov Chain Monte Carlo Stochastic Approximation Algorithm
(MCMCSAA) was developed to compute estimates of parameters with incomplete
data. In theory this algorithm guarantees convergence to the expected fixed points.
However, due to its flexibility and complexity, care needs to be taken for imple-
mentation in practice. In this paper we show that the performance of MCMCSAA
depends on many factors such as the Markov chain Monte Carlo sample size, the
step-size of the parameter update, the initial values and the choice of an approx-
imation to the Hessian matrix. Good choices of these factors are crucial to the
practical performance and our results provide practical guidelines for these choices.
We propose a new adaptive and hybrid procedure which is stable and faster while
maintaining the same theoretical properties.

7.1 Introduction

We often face incomplete data such as censored and/or truncated data in sur-
vival analysis, and unobservable latent variables in mixed effects and errors-in-
variables models. It is difficult to compute estimates of parameters such as max-
imum likelihood estimates (MLE) with incomplete data because the likelihood
usually involves intractable integrals. For example, the likelihood function of the
generalized linear mixed models (GLMM) involves an integral with respect to the
random effects which usually does not have a closed form for non-Gaussian obser-
vations. Laplace approximation may lead to large bias and inconsistent estimates
[3,7, 11]. McCulloch [13] modified the EM and the Newton—Raphson algorithms
for fitting GLMMs with integrals approximated by Monte Carlo (MC) methods. A
drawback of the MC approach is that the iterative scheme converges to a random
variable rather than to the expected fixed point. As a consequence, it is difficult
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to decide the MC sample size and a stopping rule for such an iterative procedure.
Booth and Hobert [2] pointed out these potential problems and proposed an empiri-
cal approach to increase the MC sample size. However, it is unclear if their algorithm
converges in theory.

MCMCSAA proposed by [5] (Gu and Kong, 2000, Personal communication)
for incomplete data is a major breakthrough which guarantees convergence to the
expected fixed points in theory. MCMCSAA uses stochastic approximation (SA)
to update the parameters and Markov chain Monte Carlo MCMC) to approximate
integrals at each iteration. In this article we investigate the performance of MCMC-
SAA using simulations. Our goal is to reveal some practical issues involved in the
implementation of MCMCSAA. We find that MCMCSAA can be sensitive to the
choices of (a) the initial values, (b) the MCMC sample size and step-size of the
parameter update, and (c) a matrix whose expected value approximates the Hes-
sian of the target function. We provide recommendations for implementation and
propose a hybrid algorithm which is stable and considerably faster.

We review the MCMCSAA in Sect.7.2. We conduct simulations to illustrate
potential pitfalls in the implementation of the MCMCSAA in Sect. 7.3. We propose
a hybrid algorithm in Sect. 7.4. Concluding remarks are made in Sect. 7.5.

7.2 Markov Chain Monte Carlo Stochastic Approximation
Algorithms

In this section we briefly review MCMCSAA proposed by [5] (Gu and Kong,
2000, Personal communication). See also [6, 9, 10] (Gu and Zhu, 2002, Personal
communication).

Suppose that we want to solve the following estimating equations

h(6) =0, (7.1)

where 6 is a vector of parameters and 4(6) is a vector valued function that can be

written as the expectation of a function H ((3, ¢€) with respect to a random vector &
with density function f3(é):

h(6) = E;[H(6,8)] :/H(é,é')fg(é')d?z. (7.2)

In survival analysis, for example, data might be right censored. The score function
can be written in form (7.2) [14]. Another example is a GLMM when the random
effects are considered as missing data. Integrating respect to the vector of random
effects leads to form (7.2) [5, 13, 15].

To apply the MCMCSAA, one needs to find a matrix / ((3, ¢) such that E3[1 (5 ,0)]
~ oh / 30 in the neighborhood of the solution. We consider the following three
different choices of 1 (5 ,€):
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1,(0,8) = —3H(6,¢)/06,

1(0.8) = —0H (6.8)/00 — H(0.8)H (6.)"

15(6.8) = —0H (6,8)/06 — H(6.8)H (6.8)T + Ez[H (6, 8)E;[H (6,8)]".
(7.3)

Whenever the meaning is clear we will drop the dependence on 6 and ¢ from all
I-matrices. Also for brevity we will often refer to I;, I, or I3 when we actually
mean the algorithm based on /1, I, or I3. I1 used by [1], is usually well-behaved.
For example, it is positive definite for convex target functions. Gu and Kong (2000,
Personal communication) proposed I, and claimed that it is more efficient. /3 moti-

vated by the actual derivative of h (é) with respect to 6T [12] is new. Convergence
of the algorithms are guaranteed for all three choices of I-matrices [1,5]. (Gu and
Kong, 2000, Personal communication).

Let {yx, k > 1} be a sequence of real numbers, and {my,k > 1} be a sequence
of positive integers which satisfy the following conditions:

(C1) yx <Iforallk

(€2) ZZO=1 Vi = 00

(C3) > pz,vit¢/my < oo, for some € € (0,1)
(C4) Y32 lyk/mk — Y1/ mi—1| < o0

At iteration k, a MCMC sample of size my with equilibrium distribution
f5(é) is drawn. See [4] for an overview of the Metropolis—Hastings algorithm.

Let E,(Cl), . ,E,(ka) be the MCMC sample of size my after some burn-in. The
MCMCSAA updates the parameter vector 6 and a matrix I" as follows:

e = (1= yi) D=1 + vk,

o - (7.4)
Ok = Ok—1 + I " Hy,
where
- mg )
He =Y H(Gr. )/ mr (7.5)
j=1
mpg R .
Te =Y 1018/ my. (7.6)
j=1

The I matrix in (7.6) may take any form in (7.3). I'; acts as a proxy of the Hessian
matrix and is updated as parameters. yi is the step-size of the parameter updates.
The innovation of MCMCSAA is the introduction of two sequences {yx } and {my}.

For a constant my and y; = 1, the estimate of 6 will maintain a certain amount
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of variation along iterations due to estimating H and I by averaging over ran-
dom samples. By increasing the sample size my, decreasing step-size Yk, or both,
the MCMCSAA decreases the variation in 6 as iteration increases. Gu and Kong
(2000, Personal communication) have shown that under some regularity conditions
and when my and yy satisfy conditions (C1)-(C4), ék converges to the solution of
(7.1) almost surely. The following are three combinations of my and yi that satisfy
conditions (C1)—(C4):

Gl yr = land my = mg + k?
G2 yr =1/kand mp = mg
G3 yr =1/Vkandmg =mo+k

where my is the starting MCMC sample size. We see that when y; = 1, my needs
to increase quadratically. When m, is a constant, y; needs to decrease in the order
of 1/k. G3 is a compromise between G1 and G2.

The result of (Gu and Kong, 2000, Personal communication)provides the theo-
retical justification for the MCMCSAA. However, few research has been done to
investigate its performance in practice. The simulation results in the next section
are intended to show some potential problems that may occur in practice, and to
compare different choices of parameters of the algorithm. We propose a new hybrid
procedure and conduct more simulations in Sect. 7.4.

7.3 Simulations

To implement the MCMCSAA, one needs to decide (a) the initial values éo, (b) the
MCMC sample size mj and step-size of the parameter update yg, and (c) the form
of the I-matrix. We will use simulations to show that the stability of the algorithm
and the speed of convergence depends critically on all three choices.

We use the following simple model to illustrate challenges and potential pitfalls
involved in the implementation of the MCMCSAA. Same issues arise in more com-
plex models. Consider an experiment with n = 10 binary observations from each
of m = 20 subjects. Binary observations are generate from the following simple
GLMM:

P(y; = 11b;) = exp(bi)/(1 + exp(bi)),
b N©,0), i=1,....m; j=1,....n, (7.7)

where b; are random effects. Note that there is no fixed effects in model (7.7). The
problems we are going to show in this section are usually associated with the esti-
mation of variance components such as 6, while the estimation of fixed effects is
stable and converges quickly. Thus we set them to zero to simplify the exposition.
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Let 5 = (Vi1 ¥ine YmioYmn) and b = (b1.-++ . bm). The log-
marginal distribution of  is

15(5:6) = tog [ 55616 exptlyb:6)}d, .8

where 'fil 3 (l; ; 0) is the conditional distribution of y conditional on band! 3 (l; 10) s

the log-density of b. It can be shown that under some regularity conditions,

3l (b 9)

(5, 60) (b 0) - -
y _ [ % ] _E.
_/ Sy b)db = By s =

20 20

where fEIfz is the posterior density of b given y. Thus we can apply MCMCSAA
with e = I;|)7 and H(G, I;) = dl; (l; 0)/00. It is easy to check that for model (7.7),

H(0.b) = —m/(20) + Y b?/(26%),

i=1
L(0.b) = —m/(26%) + ) _ b} /6°.
= (7.9)

i=1

m 2
1(0.b) = 1(8.b) - [—m/(w) + Zb?/(zez)} ,
I3(6,b) = 1(6, b) + (B H (6, b))%,

Since 6 is non-negative, we set 6 = 0r_ if the update 01 + v I' ,:ll:lk < 0.
We estimate El§|y*1j1(9’5) by 1:1(9,13) = ZT:I H(G,E(j)), where b are

MCMC samples. We use the single component Metropolis—Hastings algorithm to

sample from fglﬁ (l;) with the length of burn-in set to 300 [4, 8]. Specifically, at the

pth iteration of the MCMC, we update each element b? ), e b,(np ) one by one as
follows: generate X from N(bl-(p _1), 0.56;_1) as a candidate for bl-(p ) and U from
Unif(0,1); set b = X if U < min{l,7(X)/x(b" ")} and b? = pP~V
otherwise.

We simulate data with three true values of 8: 0.5, 1.0 and 2.0. Denote 6, as the
initial value and 6y r as the MLE. We compute 6,z s numerically by maximiz-
ing (7.8) using integration and optimization functions in R (www.r-project.org). We
consider the following combinations of parameters for the algorithm:

Initial values 901 0.50y1E, Ovrr and 20y £
I -matrices: I, I, and I3

yx and my: G1, G2 and G3

mo: 30 and 300
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Fig. 7.1 Four patterns of sequences divided by Oy

We have also tried to set both m and burn-in period to 10,000, the convergence
behavior remains the same. For each combination of parameters, we repeat simula-
tion 100 times. Following (Gu and Kong, 2000, Personal communication), we stop
the algorithm after 50 iterations for G1, 1,000 iterations for G2, and 250 iterations
for G3.

We first look at the convergence behavior. All iteration sequences follow one of
the following four patterns: (a) converged to the MLE, (d) approaching but not yet
converged to the MLE, (c) converges to zero, and (d) diverges to infinity. We show
a typical example for each pattern in Fig. 7.1.

The curve of sequences usually becomes smoother and smoother along the iter-
ations. However, depending on the choice of y; and mj, random variation persists
even after a large number of iterations. The following definitions allow the estimates
to have small variations around the MLE. Let 65 be average of the estimate at the
last 5 iterations. For any sequence, we define the following three states:

(S1) converged, if |(é5 — Omie)/ Opre + 1)| < 0.05

(SZ) diverged, l_f |(95 — 9MLE)/(9MLE + 1)| > 1, or |(95 — QMLE)/(QMLE + 1)| >
0.05 and |65 /6Op1e| < 0.05

(S3) not converged, otherwise

Not converged sequences (S3) consists of two classes: sequences that are converging
but are not close enough to the MLE and sequences that are diverging but are not
far enough from the MLE. Table 7.1 shows the convergence behavior. An entry i /j
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Table 7.1 Number of simulation replications that are converged or diverged

0 9() my = 30 mgy = 300
Gl G2 G3 Gl G2 G3
I 78/0 0/0 72/0 78/0 0/0 75/0
0.50m1E L 8/85 55/26 43/51 11/86 74/17 61/32
I; 38/47 53/6 70/15 50/38 80/6 75/15
I 79/0 64/0 85/0 83/0 92/0 89/0
0.5 160m1E L 5/93 55/36 32/64 21/75 84/12 62/30
I; 38/54 65/8 60/18 53/36 85/9 71/14
I 82/0 46/0 87/0 87/0 55/0 90/0
1.560m1E L 4/95 27/65 16/80 11/88 46/47 24/71
I; 34/54 63/9 50/36 50/44 83/5 71/22
I 95/0 0/0 94/0 97/0 0/0 94/0
0.50m1E I 10/85 65/22 49/44 22/77 7117 72/24
Iz 79/19 75/3 83/6 79/17 97/1 92/2
I 97/0 86/0 98/0 95/0 100/0 99/0
1 160mLE I 9/87 66/29 58/38 51/48 96/3 89/9
Iz 64/33 81/6 81/7 81/18 98/0 94/6
I 96/0 77/0 98/0 97/0 89/0 98/0
1.560m1E I 3/95 28/64 26/72 6/93 38/54 30/69
Iz 67/31 82/6 86/9 80/19 89/3 9177
I 98/0 0/0 98/0 98/0 0/0 99/0
0.50mLE L 11/88 66/24 58/40 32/66 53/42 49/49
I; 84/14 95/1 97/1 91/9 100/0 100/0
I 98/0 97/0 99/0 99/0 100/0 100/0
2 160m1E L 12/87 85/13 58/40 58/42 99/1 95/4
I; 85/15 96/0 93/5 91/8 100/0 99/1
I 98/0 87/0 99/0 100/0 98/0 100/0
1.560m1E L 8/91 35/64 19/81 5/95 16/84 12/88
I; 82/17 78/9 91/8 92/5 82/3 98/0

indicates that there are i converged sequences, j diverged sequences, and 100—i — j
not converged sequences.

It is clear that the convergence behavior depends on 6 as well as the choices of
I, yx and my. As always, a good initial value is important. /; is stable as expected:
most all sequences based on /; converged and none of them diverged. A closer look
indicates that all not converged sequences are actually converging slowly. In contrast
to /1, I and I3 are unstable with many sequences diverged, regardless of the choice
of 8y, I, yx and my. It is important to note that the problem persists even when the
initial value is set to the target value 8y . I» has more diverged sequences than /3.
Convergence also depends on the specific combination of I, I», I3 and G1, G2, G3.
For example, I, always performs best with G2. And when 6y is not close to Oy,
I, performs better with G1 than with G2.

To illustrate the cause of divergence when I, and /3 are used, for a typical
diverged case, we plot 6 and I" for the first 10 iterations in Fig.7.2. We see that



104 Y. Jiang et al.

00 — o ©
© - o)
D
Q'-_
N A\~ A\~ A\~ O A\ A\~
o —
I I I I I
2 4 6 8 10
[9)
Q'-_
o
m_
o
o~ o
L o
o O
o)
T —
I I I I I
2 4 6 8 10
iterations

Fig. 7.2 Effect of negative I" on estimate of 6. The bottom plot shows the value of I" based on /5.
The rop plot shows the estimate of € at the corresponding iteration, and the solid line represents
the MLE

from iteration 7, due to MCMC simulation error, the I" values are negative (i.e.,
the I" matrices are not positive definite). This turns 6 away from the MLE. In most
cases sequences with such problems do not recover and diverge to infinity or con-
verge to zero. Zhu and Lee [15] noticed the same problem and proposed a hybrid
algorithm.

Next, we look at the rate of convergence. In Table 7.1 we see that for all com-
binations, the algorithm based on G2 has the most sequences that are classified as
not converged. To compare G1 and G2, we show two typical sequences of estimates
based on 11 with 6y = Oy and 6y = 0.560 in Fig.7.3.

The left plot shows that /0y g are close to 1 for both G1 ad G2, which means
that both sequences settle around the MLE, but the curve based on G2 is much
smoother than the curve based on G1. The right plot shows that the sequences based
on G1 converged to the MLE in fewer than 50 iterations, whereas the sequence
based on G2 has not reached the level of the MLE after 1,000 iterations.

Plots under other settings are similar. We conclude that if the starting value is
far from the MLE, G1 reaches the level of the MLE faster than G2. On the other
hand if the estimate is already at the level of the MLE, G2 settles down much faster
than G1. These results are not surprising since for G2 the step-size of the parameter
update yy is 1/ k. Thus after only a few iteration y is very small and does not allow
a large change in 6 anymore. It takes many iterations to reach the level of the MLE.
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Fig. 7.3 Ratios between the estimates and 6, for sequences based on G1 and G2. The iteration
numbers of G1 are marked at the bottom of the x-axis and iteration numbers of G2 are marked at
the top

For G1, yi is always 1. Therefore, it can reach the level of the MLE quickly, but
remain to have random variations after the level of the MLE has been reached. G3
is a compromise between G1 and G2 (not shown).

Above simulation results illustrate advantages and disadvantages of different
combinations of the / matrices, step size yx and MCMC sample size my. These
results can be used as a guide to make an algorithm stable and efficient. Overall,
we recommend /; for stability. One may use G1 and then switch to G2 after the
sequence has reached the target level (Sect. 7.4). When I, or I3 are used, one needs
to check the properties of the estimates, for example, if the I” matrices are positive
definite. One can start with /1 and switch to I, or I3 after a number of iterations
[15]. This would combine the stability of /; in the beginning of the algorithm with
the faster convergence of I, or I3 at the end of the algorithm.

7.4 A Hybrid Algorithm

We consider /; only in this section. Based on simulations in the previous section, we
found that G1 reaches the MLE quickly and G2 reduces variation in the estimates
quickly after the sequence reached the level of the MLE. Therefore, we consider the
following class of algorithms:

Ye = k7% and mg = mo + k207 0 < <1. (7.10)

Special cases include G1 with #; = 0, G3 with #; = 0.5, and G2 with #; = 1. Note
that the sequences need to satisfy conditions (C1)—(C4).
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We would like #; to be close to zero for small k& and close to one for large k.
The idea of our procedures is to select y; and mj adaptively according to the actual
convergence behavior. There could be many adaptive schemes. We consider a simple
approach by fitting a regression line through the last K estimates of 6. If the slope
is zero, it is likely that the estimates have reached the level of the MLE and the
variation of @ around the regression line is likely due to MCMC sampling. In this
case we want to reduce the variation using a combination of y; and my similar to
G2. If the slope is not zero, i.e., the sequence is still increasing or decreasing, it
is desirable to choose yj and my such that the estimates reach the level of MLE
quickly. One such choice is G1. Let r be the correlation coefficient between the
estimate of @ at the previous K iterations and their corresponding iteration numbers,
and 7y, be the sample correlation coefficient. Zero ry corresponds to the zero slope
of the regression line. We use t-test to test hypothesis Hy : ry = 0 v.s. Hy : r # 0.

Specifically, Hy is rejected when ‘fk/‘/(l — i)/ (K — 2)‘ > to/2,K—2 Where o is

the significant level and #4/5 g—» is the 1 — «/2 percentile of the z-distribution with
K — 2 degrees of freedom. This suggests the following algorithms:

G4 Fork>K,tk=1—r]§

G5 Fork > K, tx = (1 =r)I(|fx/ /(1 =72/ (K = 2)| < ta/2,k-2)
G6  Fork > K, 1y = I(|fx/ /(1 = 72)/(K = 2)| < ta/2,k-2)

G1 is used for the first K iterations in G4, G5 and G6. To implement algorithms G4,
G5 and G6, we need to decide the number K, that is, the last K estimates to be used
to compute the correlation coefficients.

For the same simulated data we apply algorithms G4, G5 and G6 with four dif-
ferent choices of K: 10, 20, 30 and 40. We stop the hybrid algorithms after the
50th iteration which is consistent with the number used for G1 in the previous sec-
tion. Table 7.2 presents the number of converged sequences out of 100 simulation
replications. There is no diverged sequences.

Table 7.2 indicates that the convergence behavior is insensitive to the choice of
K so long it is large enough (>10). To compare the convergence rate with G1, we
plot a typical replication of hybrid algorithms with K = 20 in Fig. 7.4. We see that
the curves based on hybrid algorithms are much smoother than those based on G1.
However, it is unclear whether 50 iterations are enough for the sequences to reach
the Oy.. We need to consider stopping rules.

Different convergence criteria have been used in practice. One simple rule is
to stop the algorithm when |0x4+; — x| < 8 where § is a preassigned number.
Gu and Zhu [6] proposed a two-stage algorithm where the first stage stops at K;
where K1 = inf{K > Ko : ||Z,§:K_KO+1 %H < 81}, Ko and §; are
preassigned constants and sgn is the signum function of a real number x which
returns 1 when x > 0, 0 when x = 0 and —1 when x < 0. The second stage utilizes
the trace of 1" matrix and H matrix to decide the stopping rule. We use the variance
along iterations to measure whether the sequence is stable or not. Specifically we
consider the following two stopping rules:
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Table 7.2 Number of simulation replications that are converged

[ True value 6
0.50 1.00 2.00
G4 G5 G6 G4 G5 G6 G4 G5 G6
K =10 64 76 73 90 94 96 96 98 99
0.50y1E K =20 65 78 75 89 94 97 97 97 98
K =30 72 78 76 89 94 95 97 98 100
K =40 69 75 77 93 95 96 98 99 97
K =10 83 83 84 90 95 95 99 98 99
160y K =20 73 80 83 93 96 94 97 100 98
K =30 73 82 81 91 93 96 100 99 100
K =40 82 85 79 96 94 96 99 99 99
K =10 86 84 79 92 93 99 97 99 99
1.56y1E K =20 75 86 87 91 96 98 97 98 99
K =30 75 87 86 96 93 99 99 99 100
K =40 84 83 84 93 94 95 100 99 98
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Fig. 7.4 Ratios between the estimates and 0y ¢ for sequences based on G1, G4, G5 and G6

I At the kth iteration, stop if % < &, where 8;, 6, are predetermined

numbers and v is the sample variance of {6, ...0 }.

II Since Fk_l is an estimate of the covariance matrix of é, therefore, instead of v,
we stop if 16 —b—11
VI 8

For the same simulation settings, we apply algorithms G1-G6 using above two stop-
ping rules. For both rules, we set §; = 0.001 and 6, = 0.001. We compute k as
the average of number of iterations needed to satisfy the stopping criterion, conv. as
converged cases after 600 iterations, diff as the average of the difference between

< 82.
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Table 7.3 Convergence behavior of G1, G3, G4, G5, and G6 under stopping rule I, diff are
multiplied by 1,000 for ease of presentation

0 6o Gl G4 G5 G6 G3 |
0.5 0.50u1E k 118.59 50.12 55.02 53.53 77.40
conv. 87 68 78 78 47
diff 6.672 19.60 12.55 14.99 38.5
CPU (s) 66.12 1.34 4.38 4.67 2.64
0.5 10y k 216.40 80.38 82.44 71.86 179.42
conv. 92 82 83 84 84
diff 2.677 10.66 8.424 9.49 7.407
CPU (s) 390.82 2.71 16.49 9.80 6.77
0.5 1.5001 k 131.41 45.04 53.75 51.84 87.12
cony. 92 73 78 84 70
diff 5.881 13.90 11.46 10.16 15.23
CPU (s) 113.02 0.94 4.09 4.10 3.04
1 0.50u1E k 125.4 45.8 49.49 43.13 66
conv. 95 87 95 91 65
diff 7.588 22.13 14.15 18.86 43.98
CPU (s) 84.55 1.03 2.70 2.03 2.25
1 10y k 289.66 90.9 89.66 77.59 222.02
conv. 98 94 97 98 99
diff 3.188 12.41 9.512 11.95 10.35
CPU (s) 828.94 3.03 17.29 10.797 8.9
1 1.5001k k 156.46 90.9 56.05 49.42 88.16
cony. 98 94 95 93 89
diff 6.854 20.41 17.67 17.10 20.92
CPU (s) 199.19 1.11 4.56 3.38 3.11
2 0.50u1E k 132.87 44.82 49.33 43.01 68.16
conv. 100 97 96 98 84
diff 13.46 43.17 34.76 31.84 57.68
CPU (s) 111.41 0.93 2.92 1.88 2.31
2 10y k 272.06 96.15 90.54 79.69 239.70
conv. 99 97 100 100 94
diff 8.389 26.86 17.34 17.89 20.89
CPU (s) 720.95 3.27 14.30 11.57 9.47
2 1.501E k 162.57 49.47 55.96 47.83 80.69
conv. 100 94 97 99 94
diff 15.68 40.31 30.07 30.78 37.05
CPU (s) 195.29 1.07 4.44 2.99 2.80

estimates and the 0z among converged sequences, and CPU as the average CPU
time in seconds for one simulation replication.

Tables 7.3 and 7.4 show convergence behavior under rules I and II respectively.
We did not include the convergence behavior of G2 because G2 performs poorly
under these two stopping rules. G2 makes the algorithm stop early even though the
iteration is far away from the targeted G g.
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Table 7.4 Convergence behavior of G1, G3, G4, G5 and G6 under stopping rule II, diff are
multiplied by 1,000 for ease of presentation

0 6o Gl G4 G5 G6 G3 |
0.5 0.50u1E k 72.58 38.59 43.95 39.79 43.32
conv. 83 59 70 70 24
diff 9.562 23.15 17.69 18.51 59.47
CPU (s) 14.02 1.55 3.81 2.55 1.40
0.5 10y k 78.08 32.16 34.53 32.79 35.41
conv. 88 72 75 77 73
diff 7.743 16.12 15.23 13.35 14.72
CPU (s) 16.28 1.20 1.76 1.68 1.16
0.5 1.5001 k 68.98 32 35.08 34.32 38.89
cony. 88 74 77 76 57
diff 8.929 16.53 13.36 14.36 26.09
CPU (s) 11.97 1.18 1.85 1.77 1.27
1 0.50u1E k 71.37 33.06 38.46 36.41 38.48
conv. 96 81 91 91 41
diff 11.90 28.44 21.90 24.23 66.73
CPU (s) 13.83 1.26 2.17 1.96 1.24
1 10y k 63.86 30.2 33.91 32.68 35.98
conv. 94 87 91 89 92
diff 11.16 23.01 18.18 20.98 19.49
CPU (s) 10.07 1.12 1.68 1.61 1.17
1 1.500m1 k 65.60 30.88 31.86 33.01 36.02
cony. 98 92 91 88 84
diff 10.75 21.11 21.80 24.16 27.19
CPU (s) 12.44 1.14 1.48 1.64 1.17
2 0.50u1E k 67.01 34.21 34.19 32.87 35.21
conv. 98 94 96 97 61
diff 22.34 48.81 43.22 42.02 13.57
CPU (s) 11.25 1.28 1.66 1.62 1.14
2 10y k 65.75 29.83 29.85 27.5 32.81
conv. 100 96 96 97 95
diff 21.46 47.51 39.91 48.30 37.52
CPU (s) 10.89 1.10 1.30 1.13 1.07
2 1.501E k 68.42 29.67 32.35 30.73 33.84
conv. 100 95 99 96 99
diff 23.19 50.40 41.64 47.49 51.93
CPU (s) 12.85 1.10 1.53 1.38 1.10

First, we see that stopping rule I is much more time-consuming than stopping
rule II for G1, but not for the hybrid algorithms. And when 6y = 6Oyg, G1 with
stopping rule I is very slow to converge, the average CPU time is almost 200 times
of those for hybrid algorithms.

Overall, G1 has the highest converged cases among all the algorithms. On the
other hand, G1 is always the most time-consuming one. G3 usually has the lowest
converged cases especially when 6 is far away from the Oy g. G3 is the tradeoff



110 Y. Jiang et al.

between G1 and G2 and the sequences based on G3 is much smoother than G1. So
if we use the variation of sequences as the stopping rule, G3 may stop prematurely.
The hybrid algorithms overcome this problem by switching between G1 and G2,
so they give better converged rate than G3 even though the average CPU time for
hybrid algorithms and G3 are at the same level.

Now we look at the average difference between estimates and 6y, . Under stop-
ping rule I, when § = 0.5 and § = 1, the average difference is at 10~ level for
G1, and at 1072 level for hybrid algorithms; when 68 = 2, the average difference
is at 1072 level for both G1 and hybrid algorithms. Under stopping rule II, when
6 = 0.5, the average difference is at 1073 level for G1, and at 1072 level for hybrid
algorithms; when 6 = 1 and # = 2, the average difference is at 1072 level for both
G1 and hybrid algorithms. The average difference based on G1 are always smaller
then the hybrid algorithms. However, the CPU time for G1 is between 10 to 200
times of that for the hybrid algorithms.

7.5 Conclusions

MCMCSAA is a powerful tool to compute estimates of parameters with incomplete
data. It has attractive theoretical properties. However, due to its complexity and flex-
ibility, the implementation of MCMCSAA is far from straightforward and caution
needs to be exercised.

The MCMCSAA involves several parameters including the form of the /-matrix,
the MCMC sample size my and step-size of the parameter update y;. We have
shown that the performance of MCMCSAA depends critically on all three param-
eters. We recommend [; over I, and I3 for stability. When I, or I3 is used, one
should check the property of the H matrix to avoid divergence. There can be many
choices for the combination of the MCMC sample size my and step-size of the
parameter update yi. Different choices represent different compromise between
convergence rate and CPU time. There is no ultimate best algorithm for both pre-
cision and efficiency. Therefore, different choice should be chosen for different
purpose. In general, the proposed hybrid algorithms are stable and fast.
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Chapter 8
Stochastic Comparisons of Spacings
from Heterogeneous Samples

Subhash Kochar and Maochao Xu

Abstract In this paper we review some of the recently obtained results in the
area of stochastic comparisons of sample spacings when the observations are not
necessarily identically distributed. A few new results on necessary and sufficient
conditions for various stochastic orderings among spacings are also given. The paper
is concluded with some examples and applications.

8.1 Introduction

Spacings are of great interest in many areas of statistics, in particular, in the char-
acterizations of distributions, goodness-of-fit tests, auction theory, life testing and
reliability models. A large number of goodness-of-fit tests are based on functions of
sample spacings (see [2, 14, 15]).

Let Xy, ..., X, be n nonnegative random variables. The random variables D;., =
Xin—Xi—tpand D}, = (n—i+1)D;,i = 1,...,n, with Xo., = 0, are respec-
tively called simple spacings and normalized spacings. In the reliability context they
correspond to times elapsed between successive failures of components in a system.
In stochastic auction theory, D,., and D,., are of particular interest, which repre-
sent auction rents in buyer’s auction and reverse auction in the second-price business
auction (see [37]). It is well known that the normalized spacings of a random sample
from an exponential distribution are independent and identically distributed (i.i.d.)
random variables having the same exponential distribution. Such a characterization
may not hold for other distributions or when the observations are not independent
and identically distributed.

In many cases, the observations are independent but not identically distributed
and we call them as heterogeneous samples. The study of heterogeneous samples is
of great interest in many areas. For examples, in engineering, a complex engineering
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system is often composed of many different types of electrical components. Investi-
gating the reliability of such system is relying on heterogeneous samples, which are
the failure times of electrical components collected from experiments. Accordingly,
the topic of heterogenous samples plays an important role. However, such a study
is often challenging as the distribution theory of spacings when the observations are
not i.i.d. is often complicated. A powerful tool to investigate the stochastic proper-
ties of spacings of heterogeneous samples is stochastic orders, which is a widely
studied concept in probability and statistics.

In this paper, we review some recent results on stochastic orderings between
spacings of heterogeneous samples, and some new results and applications are
presented as well. The other interesting topic not discussed in our paper is that
of dependence among spacings. Interested readers may refer to recent papers by
[4,5,13,16,30] and references therein on this topic.

8.2 Stochastic Orders

Let X and Y be two random variables with distributions F and G, and survival
functions F = 1 — F and G = 1 — G. If X is less likely than Y to take on large
values, then, intuitively, the survival function of X will be smaller than the survival
function of Y at any fixed point. This leads to the usual stochastic order.

Definition 8.1. i( is saich be smaller than Y in the usual stochastic order, denoted
by X < Y, if F(x) < G(x), or equivalently, F(x) > G(x).

Suppose that X and Y are life lengths of two electronic components and satisfy
X <s: Y.If both components are observed to be alive at time ¢ > 0, one might con-
jecture that the residual lives would also be stochastically ordered. However, such
a result does not hold (cf. [24,26])! Hence, a stronger concept than usual stochas-
tic order is needed. The following order is motivated by the fact that the above
conjecture is false.

Definition 8.2. X is said to be smaller than Y in the hazard rate order, denoted by
X <pr Y, if

PX>x+t|X>t) <P >x+t|Y >t), forallx >0andallz.

Now, suppose that two components X and Y failed before observation time ¢ >
0.If X <y Y, is it necessarily true that the ‘inactive time’ [t — X|X < f] of X
is stochastically larger that the ‘inactive time’ [t — Y |Y < t] of Y ? The answer is
negative. The following definition is proposed to resolve this question.

Definition 8.3. X is said to be smaller than Y in the reverse hazard rate order,
denoted by X <, Y, if

Pt—X>x|X<t)>P(t—-Y >x|Y <t), forallx > 0andallzt.

An interesting order based on the mean residual life is defined as follows.
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Definition 8.4. X is said to be smaller than Y in the mean residual life order,

denoted by X <,,,; Y, if
E(X;) <E(Y;) forallt.

There are other situations, where one would like to have
[X|X € E] < [Y|Y € E]
for all possible events E. It turns out that this requirement leads to the likelihood
ratio order as defined below.
Definition 8.5. X is said to be smaller than Y in the likelihood ratio order, denoted
by X <5, Y, if
P(X >xla<X <b)<P(Y >xla<Y <b) forallxanda < b.

When the density functions of X and Y exist, X <;, Y is equivalent to

f(H)g(s) < f(s)gt) foralls <t.

The following chain of implications holds among the above univariate stochastic

orders:
Xfer=>X§th=> XfmrlY

N8 U U (8.1)
X< Y= X <Y = E(X) <E(Y).

An other order closely related to the likelihood ratio order is the joint likelihood
ordering introduced by [34].

Definition 8.6. For a bivariate random variable (X, Y), X is said to be smaller than

Y according to joint likelihood ordering, denoted by X < Y if and only if

Elp(X. V)] = E[p(Y. X)], ¢ € Gy,

where

r:{¢:¢(y’x)§¢(x’y)7 xfy}
It can be seen that
Lr:j
X1<X2 & feGy,

where f'(-,) denotes the joint density of (X1, X>).

As pointed out by [34], joint likelihood ratio ordering between the components
of a bivariate random vector may not imply likelihood ratio ordering between their
marginal distributions unless the random variables are independent, but it does
imply stochastic ordering between them, that is,

Lr:j st
X<Y=X<Y.
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A bivariate function ¢ € Gy, is called arrangement increasing (Al). Hollander
et al. [11] have studied many interesting properties of such functions, though,
apparently, they did not relate it to the notion of likelihood ratio ordering.

The above idea can be extended to compare the components of an n-dimensional

vector X = (X1,...,Xy). We define X, Zréj Zréj X, if the joint den-
sity f(x1,...,x,) of X is an arrangement increasing function. (See, [24] for the
definition of an arrangement increasing function on R”.)

In a different context, [31] studied a subclass of arrangement increasing functions
on R”, which they call as ISO* functions, as described below. Let x and y be two

vectors on R” such that ) /_,y; < >/_x; j =1,...,.n—land Y /_,yi =
> 1xi. We shall denote this partial ordering between the vectors by x < y.

Definition 8.7. A real-valued function ¢ defined on a set A C R”" is said to be
ISO* on Aif p(x) < @(y), V xKLy.

As mentioned earlier, an ISO* function is arrangement increasing but the con-
verse is not true. It is easy to see that the joint density f(x1,x2) of a bivariate
random vector (X1, X3) is ISO* if and only if the conditional density of X, given
X1 + X, =t is monotonically increasing for each fixed .

The usual likelihood ratio order has the following multivariate version.

Definition 8.8. Let X = (X1, X5,...,X,) and Y = (Y1,Y2,...,Y,) be two n-
dimensional random vectors with absolutely continuous [or discrete] distribution
functions and let f and g denote their continuous [or discrete] density functions,
respectively. Suppose that

S®gy) = fxAy)gxVy)

for every x and y. Then X is said to be smaller than Y in the multivariate likelihood
ratio order (denoted as X <;, Y).

It is known that multivariate likelihood ratio order implies component-wise like-
lihood ratio order. For more details on the univariate and multivariate stochastic
orders (see [26,33]).

A basic concept for comparing variability among distributions is that of disper-
sive ordering as defined below.

Definition 8.9. X is said to be less dispersed than Y, denoted by X <, Y, if
F'B) -F ') =G '(B) -G ()

forall0 < @ < B < 1, where F~! and G~! denote their corresponding right
continuous inverses.

For more discussion, one may refer to Sect. 3.B of [33].



8 Stochastic Comparisons of Spacings 117

A weaker order called the right spread order in [9], or excess wealth order in [32]
has also been proposed to compare the variabilities of two distributions.

Definition 8.10. X is said to be less right spread than Y, denoted by X <ggs Y, if

o0 o o0 o
/ F(x)dx < / G(x)dx, forall0<p <I1.
F~1(p) G~ l(p)

It is known that

X <aip Y = X <ps Y = Var(X) < Var(Y).

8.3 Spacings

As mentioned earlier, the normalized spacings of a random sample from an expo-
nential distribution are i.i.d. random variables having the same exponential distribu-
tion. Many authors have studied the stochastic properties of spacings from restricted
families of distributions. Barlow and Proschan [3] proved that if X;,..., X, isa
random sample from a decreasing failure rate (DFR) distribution, then the succes-
sive normalized spacings are stochastically increasing. Kochar and Kirmani [20]
strengthened this result from stochastic ordering to hazard rate ordering, that is, for
i=1,....,.n—1,

Dl*n Zhr D;k+1:n' (82)

The corresponding problem when the random variables are not identically dis-
tributed has also been studied by many researchers. For a review of this topic see
[19]. In the following, we review some important results obtained recently on this
topic and also give some new results.

First, let us introduce the proportional hazard rates (PHR) model, which will be
discussed extensively in the paper. Independent random variables X1, X2, ..., X»
are said to follow PHR model if fori = 1,2, ..., n, the survival function of X; can
be expressed as,

Fi(x) = [F()), ford; >0, (8.3)

where F(x) is the survival function of some random variable X . If r(¢) denotes the
hazard rate corresponding to the baseline distribution F, then the hazard rate of X;
isA;r(t),i =1,2,...,n. We can equivalently express (8.3) as

Fi(x) = e MR™ i — 12 . .n
where R(x) = f_xoo r(t)dt, is the cumulative hazard rate of X. The PHR model

includes many well known distributions, such as exponential, Weibull, Pareto and
Lomax, etc.
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8.3.1 One-Sample Problem

Pledger and Proschan [29] considered the problem of stochastically comparing the
spacings of heterogeneous samples in the PHR model.

Theorem 8.1. ile, ..., Xp are independent random variables with X; having sur-
vival function F(t) = e RO i = 1,... n, where R(t) is a concave (i.e., F is
DFR) and differentiable hazard function, then

DY, <ot D}, for i<,

Kochar and Kirmani [20] proved that the joint density of the normalized spacings
is ISO* when the joint density of the parent observations is convex. This will hold,
in particular, when the X;’s are independent (but not necessarily identical) with
log-convex densities.

Theorem 8.2. Let the joint density fx(x1,...,xn) of X = (X1,..., Xy) be convex.
Then the joint density of D = (D1, ..., Dpy) is ISO*.

This result immediately leads to the following interesting result.

Theorem 8.3. Let X1, X»,...,Xn be independent random variables with log-
convex densities. Then

" Lr:.j " Lr.j Lr.j "
Dl:n = DZ:n = ... = Dn:n'

If a density is log-convex, it is DFR, but the converse is not true. Hence, Theo-
rem 8.3 establishes a stronger ordering between the normalized spacings than does
Theorem 8.1 under a stronger condition on the parent distributions. A related paper
on this topic is by [27].

One may wonder whether under the condition of Theorem 8.3 the result can be
extended from joint likelihood ratio order to likelihood ratio order? The answer is
negative as illustrated by a counterexample in [25].

Let Xy,..., X, be independent exponential random variables with X; having
failure rate A; fori = 1,...,n. Hu et al. [12] proved the following result.

Theorem 8.4. If A, +1 < [>]Ar fork =1,...,n, then
Dy =i Dpyinst

and
Dl:n <Ir D2:n+1[D2:n+1 <Ir D2:n]-

IfA;i + Aj > Ay for all distinct i, j and k then

Dn—l:n <Ir Dn:n, Dn:n+1 <Ir Dn:n~
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They also showed that D,.3 <;, D3.3 forall A;’s, and
Dy <ty Doy, n>2.

Observing (8.2), [21] posed the following interesting question. Let X1, ..., X,
be independent exponential random variables with possibly different scale parame-
ters. Will it hold that

D}, <pr Dy, i=1...n-1? (8.4)
They proved their conjecture for n = 3, but the general case still remains an open
problem. This topic has been extensively studied by [6, 18, 35, 36] in single-outlier
or multiple-outlier exponential models.

8.3.2 Two-Samples Problem

Pledger and Proschan [29] considered the problem of stochastically comparing
the spacings of nonidentical independent exponential random variables with those
corresponding to stochastically comparable independent and identically distributed
exponential random variables.

Theorem 8.5. If X1,..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n,andYy,..., Y, isa mn_dom sample of size
n from an exponential distribution with common hazard rate AL = Y '_, Ai/n,
then

H*n Sst Dl*n

1

where H, = (n —i + 1)(Yj;n — Yi—1:) for i = 1,...,n are the normalized
spacings of Y;’s, and Yo., = 0.

Kochar and Korwar [21] strengthened and extended this result from stochastic
ordering to likelihood ratio ordering and dispersive ordering.

Theorem 8.6. If X1,..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n,andYy,..., Y, isa mn_dom sample of size
n from an exponential distribution with common hazard rate AL = Y '_, Ai/n,
then

H =ir Dy,

* *
in in and Hi:n Sdisp D;

e =100

Kochar and Rojo [22] further strengthened Theorem 8.6 to multivariate likeli-
hood ratio order.

Theorem 8.7. If X,,..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n,and Y1,...,Y, isa ran_dom sample of size
n from an exponential distribution with common hazard rate AL = Y i_, Ai/n,
then,
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(Hln""’HrT:n) =ir (Dik:n“"’D;::n)

As a consequence, [22] pointed out that
Yim —Yin <st Xjin — Xizp forall 1=<i<j=<n.

Let Xy,..., X, be independent exponential random variables with X; having
hazardrate A;,i = 1,...,n,and Y7, ..., Y, be a random sample of size n from an
exponential distribution with common hazard rate A. One natural question is to find
sufficient and necessary conditions for comparing simple sample spacings of Dy,
from X;’s and Hg., from Y;’s. Kochar and Korwar [21] proved that the distribution
function of D, fori = 2,...,n is a mixture of independent exponential random
variables Wlth the density functlon

[Ti=y A (Z;lfk Ar ) exp { - Z’Jl'=k Ars

)

* X) =
ka:n() gﬂ?zlz'}:ikrj n—i+1 n—i-+1

where r extends over all of the permutations of {1, 2, ..., n}. Hence, the distribution
of Dy, could be represented as

FDk:n (X) = Z ij)L:j (X),

JEr
where j denotes a permutation of (A1, ..., A,) belonging to r and

P = [T7=y A
SR EEDY R

and

* Z;l'=k Ar/

K™=k + 12
and F’ Ay, means an exponential distribution with hazard rate )&*
Note that the distribution function of Hg., is

Ax
FHy, () = Faj—k+1)(x) = 1 — eXp{_n —k+ 1%'

The following results provide sufficient and necessary conditions for stochasti-
cally comparing Dg., and Hg., according to likelihood ratio and reverse hazard rate
orderings.

Theorem 8.8. Let X1, ..., X, be independent exponential random variables with
X; having hazardrate A;, i = 1,...,n, and Y1, ..., Y, be a random sample of size
n from an exponential distribution with common hazard rate A. Then, for k > 2,

(a) Hi:n <1y Dg:p or
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(b) Hk:n =rh Dk:n
if and only if

H?= A,‘ n 2
1 2 m (Zj=k Arj)

>
Tn—k+1 vy Mgk oy
> H?:IZTEI-:,- i, D=k Ar;

Proof. The necessary and sufficient condition for likelihood ratio order between
Hpy., and Dy, follows from Lemma 3.1 of [40]. It remains to prove that the
condition on A is a necessary condition for the reverse hazard rate order.

Note that the reverse hazard rate of Dy, for k > 2, by Taylor’s expansion around
zero, is

P (x) = Ljer Piiy exp{_kl}ijx}
) ZjErpj(l —CXP{—AkJ-X})
_ Zjer pjlltj(l - A]tjx +0(x))
Zjerpj/x]tjx +o(x)
_ Zjer pjkltj - Zjer pj(/lzj')zx + o(x)
Zjapj/\zjx + o(x)

Similarly, fori > 2,

A—=A%x/(n—k 4+ 1)+ o(x)

PHin () = Ax + o(x)

Then
FHy., (X) < Fpy., (X)
implies
2
Zjer pj (M:j)
A>(m—k+1)————7—.
Zjer pJAkj

The result follows immediately. O

Analogously, we have an equivalent necessary and sufficient condition for hazard
rate order, dispersive order and stochastic order between Dy, and Hy.,,.

Theorem 8.9. Let X1, ..., X, be independent exponential random variables with
X; having hazard rate A;, i = 1,...,n,and Y1, ...,Y, be a random sample of size
n from an exponential distribution with common hazard rate A. Then, for k > 2,

(a) Hi:n <pr Di:n or
(b) Hi:n =disp Dy, or
(¢c) Hi:p <st Di:n
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if and only if

i=1
zn_k+1zn—1211 Zkr/

rjjk

Proof. Using Lemma 2.1 in [28], it is easy to show the condition on A is a neces-
sary and sufficient condition for hazard rate order between Dy., and Hy.,. Since
Hy.,, is an exponential random variable, according to Theorem 3.B.20 of [33], (a)
implies (b). By Theorem 3.B.13 there, (b) implies (c). Hence, it is enough to show
the condition on A is the necessary condition for the stochastic order. Using Taylor’s
expansion around zero for the distribution function, for k > 2,

Fpi,,(X) =Y pi(Af;x + 0(x)),

JEr
and
FHk:n (X) = mx + O(X).
So,
FHk:n (X) 2 FDk:n (X)
implies,
A=m—k+1)Y piAr.
jer

Hence, the required result follows. O

Below is another result in the same direction.

Theorem 8.10. Let X1, ..., X, be independent exponential random variables with
X; having hazard rate A;, i = 1,...,n, and Y1, ..., Y, be a random sample of size
n from an exponential distribution with common hazard rate A. Then, for k > 2,

(a) Hi:n <mri D or
(b) Hx.y <Rrs Dp:p or
(¢) EHy.p < EDpg.y

if and only if
1 1
Sk T
n— i=1
Z Z/ k)W H[ IZ/ IA”‘

Proof. From Lemma 2.6 of [38], it is seen that the condition on A is necessary and
sufficient for the mean residual life order. By Theorem 3.C.6 of [33], (a) implies (b).
It is easy to see that (b) implies (c). Hence, it is enough to show the condition on A
is a necessary condition for (c). Note that

E(Dk:n) Z A* ’
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and
n—k+1

1 )

the result follows immediately. O

E(Hk:n) =

Example 8.1. Suppose X1, X», X3 are independent exponential random variables
with parameters A1, A5, A3, respectively; and Y7, Y5, Y3 are i.i.d. exponential ran-
dom variables with parameter A. Then, from Theorem 8.9,

Y33 — Y2:3 <pr X33 — X23

if and only if

L >, o ikl 1 1t 1
= ot A Ut As At As M+ )

From Theorem 8.8,
Y33 — Ya:3 <jp X3:3 — X333

if and only if

A>A_3( LR SR )“
- lr_z Ar + A3 A1+ A3 AL+ A '

From Theorem 8.10
Y3:3 — Y2:3 <gps X33 — Xa:3

if and only if

AzARs=[ A1Aaks ( A2+ A2 A2 4 A2

A2z 4243 N2 +43)A325 (A +A3)A7A5
A2+ 22 )r
2T ).
(A2 + A)A3A3
It is worth noting that from (8.1), it follows that
Alr = Anr = ARS-

For example, let
A =1, =213 =3.

Then,
Ay = 1.91489 > Ap, = 1.56667 > Agps = 1.30435.
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8.4 Sample Range

Sample range is one of the criteria for comparing variabilities among distributions
and hence it is important to study its stochastic properties.
Kochar and Rojo [22] proved the following result.

Theorem 8.11. If X1, ..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n,and Y1,..., Y, is aﬁandom sample of size
n from an exponential distribution with common hazard rate . =y ¢ _; A; /n, then,

Yn:n - Yl:n Sst Xn:n - Xl:n-
The next theorem due to [17] improves upon this result.

Theorem 8.12. If X1, ..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n, and Y1, ..., Yy is a random sample of size
n from an exponential distribution with common hazard rate A = (I, )k,-)l/n,
then,

Yn:n - Yl:n Sst Xn:n - Xl:n-

Recently, [39] obtained a necessary and sufficient condition for stochastically
comparing sample ranges from heterogeneous and homogeneous exponential
samples.

Theorem 8.13. If X1, ..., X, are independent exponential random variables with
X; having hazard rate A;, i = 1,...,n, and Yy, ...,Yy is a random sample of size
n from an exponential distribution with common hazard rate A, then,

N l—[ﬂ A 1/(n—1)
Yn:n_Yl:n Sst Xn:n_Xl:n@/\ZAZ(%) ’

where X = Y1_, Ai/n.

Note that A < 5&, which improves upon the results of Theorem 8.12.
Kochar and Xu [23] partially strengthened Theorem 8.11 from stochastic order
to reverse hazard rate order.

Theorem 8.14. Let X1, ..., X, be independent exponential random variables with
X; having hazard rate A;, i = 1,...,n. Let Yy1,...,Y, be alandom sample of size
n from an exponential distribution with common hazard rate . =Y, A; /n. Then

Yn:n - Yl:n =rh Xn:n - Xl:n-

They also proved the following theorem for the PHR model.
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Theorem 8.15. Let X1,..., X, be independent random variables with X; having
—=Ai . .
survival function F°, i = 1,...,n. Letin, ..., Yy be a random sample with

. . e . =A -
common population survival distribution F~, where A = Z;’:l Ai/n, then
Yn:n - Yl:n st Xn:n - Xl:n-

More recently, [10] proved the following result.

Theorem 8.16. Let X1, ..., X, be independent exponential random variables with
X; having hazard rate A;j, i = 1,...,n, and Y1,...,Y, be a random sample of
size n from an exponential distribution with common hazard rate . = 3 71—, A; /n.
Let ¥y (x) and 7y (x) be reversed hazard rates of Xp.y — X1:n and Y.y — Y1,
respectively. Then

Fx (x)

7y (x)

is increasing.

The following results follow as consequences as shown in that paper.

Corollary 8.1. Let X1,..., X, be independent exponential random variables with
X; having hazard rate A;, i = 1,...,n. LetYy,...,Y, bea }Emdom sample of size
n from an exponential distribution with common hazard rate A, then,

Yn:n - Yl:n =Ir Xn:n - Xl:m

and
Yn:n - Yl:n fdisp Xn:n - Xl:n-

8.5 Applications

8.5.1 Type-I1I Censoring

If n items are put on life test and the test terminated at the time of rth failure, then

r r
— 7 — *
- 2 : - RO —I - E : in
Irn (n 1+ 1)(X,n Xl' 1,,) D

i=1 i=1

represents the total time on test (TTT), which is a well-known concept in statistical
reliability. As pointed out in [8], if those component lifetimes are i.i.d. exponential
with mean 6 or equivalently the failure rate A = 1/6, the best estimator (UMVUE)
of 0 is
h

pt

0 =

It is well-known that (cf., [8]),
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2Trn L,
— X2r>

r

where 3, denotes a chi-square distribution with 2r degrees of freedom. Based on
this result, many estimation and testing statistical procedures have been established.
However, if the components are actually from different exponential distributions
with failure rates )LT, ..., Ay, then it follows from Theorem 8.7 and Theorem 6.E.8
of [33] that ) )

0% >4 6,

where . .
é* — Zi=1 D itn
r
and A = Z?:l )L?‘ /n. This means that based on TTT statistic, one would overesti-
mate the average lifetime of components using heterogeneous exponential compo-
nents.

8.5.2 Reliability

An n-component system that fails if and only if at least k of the n components fail is
called a k-out-of-n: F system. The lifetime of such a system could be represented as
Xi:n- The k-out-of-n system structure is a very popular type of redundancy in fault
tolerant systems with wide applications in industrial and military systems. For two
different systems say a k-out-of-n: F system and a k + 1-out-of-n: F system, the
engineer may be interested in the additional lifetime Xy 1., — X, for the system
design and the cost purpose. Due to the complicated distribution form, one may
provide a sharp bound on the survival function of this based on Theorem 8.9.

For example, a plane has four engines, and a minimum of three engines are
required for the plane work. Hence, this plane is a 2-out-of-4: F system. If the
engineer is planning to improve the system to the 3-out-of-4: F system, he/she has
to consider the cost and the reliability of improvement simultaneously. Hence, it
is important to estimate the survival probability and the mean additional lifetime
X3:4 — X2:4. Theorem 8.9 provides a lower bound for the survival function of the
additional lifetime. Let (A1, A2, A3, A4) = (1,2, 2.5, 3), then,

P(X3:4 — Xo:4 > x) > e 097%,
Using Theorem 8.10, the following sharp lower bound could be established,
E(X3:4 — X5:4) > 1.0879.

In the following table, we list the lower bounds on A’s for various partial orders
to hold in Theorems 8.8-8.10.
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A Ir/th hr/ disp/st mrl/RS/E
Dj.4 2.060897 2.039216 2.019271
D34 1988454 1909626 1.838388
D4y 2.009722  1.674501  1.382051

The values above are comparable to the arithmetic mean 2.125, geometric mean
1.96799 and harmonic mean 1.791045 of A;’s. It is interesting to note that all of
the values are less than the arithmetic mean, which coincides with the condition in
Theorem 8.7.

8.5.3 Dependence Orderings Among Order Statistics

Recently [1] proved that, in a very general sense, the dependence between pairs of
order statistics decreases as the indices of the order statistics draw apart when the
observations are independent and identically distributed. Their proofs make use of
the various results on stochastic orderings between spacings and order statistics in a
nice way.

Let Xi,..., X, be independent continuous random variables with X; having

survival function fxi, i =1,...,n,and Yq,...,Y, be ii.d. continuous random
variables. Dolati et al. [7] used the concept of more RTI (right-tail increasing)
dependence order to investigate the relative dependence between the extreme order
statistics. They showed that

(Xn:n|X1:n) <r11 Ynen|Y1on).

They wondered whether the result can be extended from more RTI dependence
order to more SI (stochastic increasing) dependence order which is a stronger depen-
dence order. Genest et al. [10] have successively made use of Corollary 8.1 to solve
this problem by proving

(Xnn|X1:n) <st Ynn|Y1:n).
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Chapter 9
The Distributions of the Peak to Average and
Peak to Sum Ratios Under Exponentiality

Tomasz J. Kozubowski, Anna K. Panorska, and Fares Qeadan

Abstract Let Eq, E,, ..., Exn be independent and identically distributed exponen-
tial random variables, and let Y = \/lN:1 E;iand S = Z;N=1 E; be their maximum
and sum, respectively. We review distributional properties of the peak to sum and
peak to average ratios, R = Y/S and R = Y/(S/N), respectively, with determin-
istic N, and provide an extension to the case where N is itself a random variable,
independent of the { £;}. Our results include explicit formulas for the relevant den-
sity and distribution functions, which apply to any distribution of N, as well as a
particular example with geometrically distributed N. An example from climatology
shows modeling potential of these models.

9.1 Introduction

Let £, E», ... be independent and identically distributed (IID) exponential random
variables with the probability density function (PDF)

f(x) = BeP*, x>0, 9.1)

and let N be a discrete random variable taking values in the set N = {1,2,3,...}
of positive integers, independent of the {E;}. Let X = ZlNzl E; be the sum and
Y= \/fv=1 E; be the maximum (peak) of the random number N of the { E;}. In this
paper we study distributions of the following two ratios: the peak to sum ratio

Y N E
R=_ — \/11;—1 9.2)
X Yi=1 Ei
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and the peak to average ratio

N
Y _ \/i=1Ei

R= = )
N
X/N %Zi:lEi

(9.3)

While our results are valid for any distribution of N, we concentrate on the particular
case where N is a geometric random variable N ~ Geo(p) with the PDF

m=P(N=n)=pl-p"', neN (9.4)

Let us note that when the number of the {E£;} is a random variable, there is no
simple relation between the distributions of the two ratios R and Ié, which is in
contrast with the case when N is deterministic.

The distributions of the rations have been studied for many years, because of
their importance in various applications, especially in engineering, communications,
computer engineering, scheduling, and insurance (see, e.g., [1,6, 17,21] and the
references therein). Perhaps the most important applications of R and R are in engi-
neering, where peak to average power ratio typically needs to be minimized, see
[21] and the references therein. However, very little is known about the exact distri-
butions of the ratios with general distribution of the { £ ; } and either deterministic or
random N, except for the case of exponential distribution and deterministic N, for
which explicit formulas are available for the PDF as well as the CDF. These results,
which actually go back to [8], appeared in the context of testing for outliers in expo-
nential samples (see [12, 15]), where the ratios of order statistics to their sum are
in common use. However, these results are not widely known. In one of the earliest
works, the authors [17] derive the exact distribution of the peak to median ratio,
arguing that using median rather than mean allows for standard derivation as both
the peak (maximum) and the median are order statistics. There are also numerous
works dealing with the asymptotic results for the ratios, as well as for the distribu-
tion of the vector (X, Y), none of which present the explicit exponential case (see,
e.g.,[2,4-6,9,10,16,18]). We hope that our brief summary of the deterministic case
with exponentially distributed { E; } will be helpful in this regard.

We consider the ratios (9.2) and (9.3) with both deterministic and random N.
We shall denote the distribution of R by PTSRn or PTSRN (peak to sum ratio),
according to whether the number of terms is deterministic or random. Similarly, we
shall use the notation PTARn or PTARN (peak to average ratio) for the distribution
of R. We first review the exponential case with deterministic number of terms in
Sect. 9.2. In Sect. 9.3 we present new general results for random N. A special case
with geometric N is treated in Sect. 9.4, while a practical illustrative data example
from climatology appears in Sect.9.5. Technical derivations are deferred to the last
section.
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9.2 Models with Deterministic Number of Terms

Here we review basic results related to the ratios R and R with deterministic number
of terms, including PDF and CDF for both ratios.

9.2.1 Peak to Sum Ratio

Let R have a PTSRn distribution defined by (9.2), where N = n is non-random
and Eq, E,, ... E, are IID with exponential distribution (9.1). Clearly, whenn = 1,
the distribution of R is a point mass at 1 with probability one. One way to obtain
the PDF of R = Y /X for general n > 2, is via the joint distribution of the sum
X = Z:’-zl E; and the maximum ¥ = \/lN:1 E; of 1ID exponentials, recently
derived in [20]. A standard transformation R = Y /X, S = X leads to the joint
PDF of (R, S), which actually turns out to be the product of the marginal PDFs of
R and S, showing the independence of these quantities — which is a well-known
property of exponential samples — as well as the exact form of the PDF of R, shown
below. The support of the distribution of R is the interval [1/n, 1], and for each
t € [1/n,1], the PDF of R is

YK a1 =512 A <r<i k=12,...n-1

ha(1) = k¥l = ©.5)
otherwise,
where
n—1 s+1
ay(s) =nn-—1) o1 (=D, (9.6)
Subsequent integration of the PDF produces the CDF, which turns out to be
0, t<1/n,
Hy(t) = {55 ()10 =—so 1, Ao <e<bik=12,...n-1,
1 t>1.
9.7)

Remark 9.1. Ttis worth noting that the quantity ay, (s) in (9.2) arises as the likelihood
ratio test statistic for an upper outlier in exponential samples (see, e.g., [7, 11-13,
15]), and its distribution was derived in this context in [15]. This was accomplished
by first establishing the recursive relation

hn(0) = nfipr(6) Hy (1#_1) Crelin 1), 9:8)
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between the PDF and the CDF of R, where

I(a+b)

a—1 b—1
F(a)F(b)t 1-0°""", telo,1], 9.9)

Jap(t) =

is the PDF of a beta distribution with parameters a, b € (0, 1). Repeated application
of (9.8) leads to the formula (9.7), which was actually derived earlier in [8] via a
rather heuristic arguments. Differentiation of (9.7) produces the PDF (9.5). Further,
as noted in [15], similar considerations apply to the ratio connected with random
samples from a gamma distribution with shape parameter o > 0, in which case the
recursive relation (9.8) still holds with f7 ,_ replaced by fy o(n—1)-

Remark 9.2. Notice that the tail probability, P(R > t), takes a particularly simple
form for ¢ € (1/2, 1), since we have P(R > t) = n(1 — )"~ 1.

Remark 9.3. Observe that when n = 2, the ratio R has a uniform distribution on
its supporting interval [1/2, 1], while for n = 3 we get a triangular distribution on
the interval [1/3, 1] with a unique mode m3 = 1/2 (see Fig.9.1). It can be shown
by elementary calculations that for n > 4 the densities (9.5) are differentiable on
the interval (1/n, 1), with the values of zero at the endpoints. It also appears that all
densities are unimodal with the unique mode in the interval [1/n, 1/2], which can
be proven by straightforward algebra for the cases n = 4, and 5, where the modes
are mg = 5/11 and ms = (30 — +/32)/62, respectively.

0 1 I
0 1/8/71/6 1/5  1/4 1/3 1/2 1
r

Fig. 9.1 The densities (9.5) of the ratio (9.2) forn = 2,3,..., 8
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9.2.2 Peak to Average Ratio

The CDF and PDF of the peak to average ratio R defined in (9.3) with N = n

(denoted by PTARn) are easily derived from those of PTSRn via the relation R 4
nR. The PDF of R is given by g, (t) = (1/n)h,(t/n), where h,(x) is the PDF of
R defined in (9.5). Note, that this PDF is nonzero for ¢+ > 1. In turn, the CDF of
PTARn is G, (¢t) = H,(t/n), where H,(x) is the CDF of PTSRn given above.

9.3 Models with Random Number of Terms

In this section, we consider a general case, were N is an integer-valued random
variable supported on the set N, with

P(N =n)=p,, neN, (9.10)

while in the following section we present a particular example where N has a
geometric distribution.

9.3.1 Peak to Sum Ratio

To obtain the CDF of the PTSRN distribution, we use a standard conditioning argu-
ment. First, observe that the probability P(R < x) is zero whenever x < 0 and one
if x > 1. Second, for any 0 < x < 1, we have

P(R<x)=) P(R=x|N=nP(N =n) =) Hny(x)pn,
n=1

n=1

with H, and p, given by (9.7) and (9.10), respectively. Assuming that 1/(k + 1) <
x < 1/k with some k € N and taking into account (9.7), after some algebra we
obtain

k %)
P(R<x)=)» (-1)° Y (Z)(l —sx)" 'pp +P(N >k +1). (9.11)
s=1 n=k+1

Note that the function (9.11) is continuous in x on the interval (0, 1), with the one-
sided limits of

x—>0"

lim P(R <x) =0, lim+ P(R<x)=1-p;.
x—>1
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The CDF of R is continuous on R except at x = 1, where there is a jump of p;.
Thus, as expected, the distribution of peak to sum ratio R is a mixture of a point
mass at 1 (with probability p;) and a continuous distribution on the interval (0, 1)
(with probability 1 — p;), with the latter given by the CDF F'(¢):

0, t <1,
F(ty={F*(1), ;47 <t<¢. keN (9.12)
1, t>1,
where
1 k > n
F*@) = 1 Z(—l)s Z ( )(l—st)"_lpn+]P’(N >k+1)
N s=1 n=k+1 §

By taking the derivative, we obtain the density of the continuous part,

k 0
_ 1 _1)\s+1 n _ _ n—
f) = =7 s§=1S( 1)*+ n=§k+1 (S)(n Ds(1=st)"2py. (9.13)

for kl? <1 < ¢,k € N. The following result summarizes this discussion.

Theorem 9.1. The CDF of R defined by (9.2) is given by
G@t)=(0—=p)F@)+ pilie(?), t €R, (9.14)
with F given by (9.12), so that
RLT+0-Dx, (9.15)

where X has the CDF (9.12) and I is an indicator variable with P(I = 1) = py,
independent of X .

9.3.2 Peak to Average Ratio

We now turn to a derivation of the PTARN distribution of (9.3). Proceeding again
with a standard conditioning argument, we obtain

P(R<x)=) P(R<xIN=mPW\N =n) =} Hu(x/n)pn.

n=1 n=1
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with H, and p, given by (9.7) and (9.10), respectively. Since H,(u) is zero for
u < 1/n, the probability ]P’(R < x) is zero whenever x < 1. Assume that x > 1,
with [x] =m € N, sothatm < x < m + 1. Since H,(u) = 1 whenever u > 1, we
have H,(x/n) = 1 whenever n < m = [x]. On the other hand, for n > m, we have
1/(k +1) <x/n <1/k with k = [n/x], so that in view of (9.7) we obtain

[n/x]
Ha(x/m) = Y (Z)(—nm —sx/ny' 7t 41

s=1

This leads to the following expression for the CDF of R:

0, 1,

G(x) = oy *=b 9.16)
Zn [x]+1 Pn ( )( DS(1—sx/n)" ' x=>1.

Note that G(7) is continuous except for a jump of p; at x = 1, since R = 1

whenever N = 1, which occurs with probability p;. Thus, the distribution of Risa
mixture of a point mass at 1 and a continuous distribution on (1, 00), the latter given
by the CDF

F(x) 0 x <1,

X) =
lpl{l_p1+z x]+1pn2n/x]()( D*(1 —sx/n)"~ 1} x> 1.
9.17)

We summarize this discussion in the following result.

Theorem 9.2. The CDF ofﬁ defined by (9.3) is given by
G(t)=(0—=p)F(@)+ pilie(?), t €R, (9.18)
with F given by (9.17), so that
RL7+(1-Dx, (9.19)

where X has the CDF (9.17) and I is an indicator variable with P(I = 1) = py,
independent of X .

9.4 Geometric Example

In this section, we illustrate our general results with an example of geometrically
distributed N with PDF given by (9.4). Let us start with the PTSRN distribution,
denoted by PTSRN(p). By replacing the quantity p, in (9.12) with the geometric
PDF (9.4), we obtain the following expression for the CDF of the random variable
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X of Theorem 9.1:

k 00
ny . a1 k-1 1 l
F(t)— g n%—l(s)(l Q)"+ (1=p), k+1§t<k,keN,
(9.20)
where
q=gqs(t;p)=1—(10—-p)(1—1s) € (0,1). 9.21)

The following result provides an explicit expression for the infinite sum in (9.20).

Lemma 9.1. Foranyq € (0,1),k € N, and s € {1,2,...,k} we have
) — k+1
1—¢q)*! —1
> (acar =2 (" ag 02
s g5t s
n=k+1 n=s+1

This leads to the result below.

Theorem 9.3. Let R ~ PTSRN(p). Then the CDF of R is given by
Gt)=(0—=p)F(t) + plo)(), t €R, (9.23)

where F(1) = 0fort <0, F(1) = 1fort = 1, while for 55 <t < .k €N,

k q)s 1 k+1 n—1
F)=— Z 1>S[ s -y ( s )(1—q>"—2}+(1—p)"—1,

n=s+1
(9.24)

with g = qs(x; p) given by (9.21).

Remark 9.4. As before, the random variable R admits the mixture representation
(9.15), where the continuous variable X has the CDF F above. Moreover, by taking
the derivative, we find the PDF of X to be

[(s +1-2¢9)(1—¢q)"2 ©9.25)

k
f)y=p Zs(—l)s+1 o

k+1
- ( )(n—le— )~ 3}

n=s+1

for kl? <t< %, k € N, and zero otherwise.

Finally, we consider the peak-to-average ratio (9.3) with geometrically dis-
tributed N, which distribution shall be denoted by PTARN(p). When we replace
the quantity p, in (9.17) with the geometric PDF (9.4), we obtain the following
expression for the CDF of the random variable X of Theorem 9.2:
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Flx) = 0, x <1,
L4 125 S g Se () D1 = p (1 —sx/m ! x : i'@

In this case the mixture representation (9.19) still holds with P(/ = 1) = p and X
having a continuous distribution on (1, co) given by the CDF (9.26).

9.5 An Illustrative Data Example

We illustrate the possible applications of the models for the ratio of the maximum
to the total of IID exponentials with an example from climatology. Understanding
the distribution of the ratio of maximum to the total precipitation over a time period
helps with water management and flood control (see e.g., [19,22,23], or [24] and
the references therein). The issue of flooding is especially important to study in the
“low countries”, such as the Netherlands. The latter is a country with about 25%
of its area below the mean sea level, making 56% of its population living on the
floodplains (see [25]) and 70% of the country gross domestic product produced in
these low areas. Through the Netherlands’ history, countless people have lost their
lives and homes to floods from the North Sea or the rivers that could not be held by
the flood defenses. The most recent flood events in the Netherlands were the 1993
and 1995 floods and the disastrous North Sea Flood of 1953 when 1,836 people
were killed (see [3]).

For this illustration, we chose precipitation data from two stations nearby a mas-
sive river delta in the southwestern Netherlands, west from Antwerp: station 745
in Axel and station 770 in Westdorpe. Axel has data records from 1906-1995 and
Westdorpe from 1995-2008. The two towns are about 4 miles apart. The first station
is located at 51°17’ latitude, 3°55’ longitude, and 2.1 (m) elevation, while the West-
dorpe station is located at 51°13’ latitude, 3°51’ longitude, and 1.5 (m) elevation.
The precipitation is measured at 08.00 UT, as a 24 h total (from 08.00 to 08.00 UT),
see "http://www.knmi.nl/"for more details. The precipitation measurement
unit is 0.1 mm. For modeling, the data consisted of 4,197 weekly precipitation totals
(from 01-01-1928 to 06-07-2008). Each four consecutive weeks were treated as
1 month. One month with zero total precipitation was excluded from analysis. We
had 1,049 months in the data set . For each month two variables were computed:
monthly total precipitation and monthly maximum precipitation. Monthly total was
the sum of the four weekly totals and monthly maximum was the largest weekly
total for the 4 weeks in the month. The data were then converted to the ratios of the
maximum to the total, resulting in 1,049 values.

We fit the distribution PTSRn with n = 4 to the precipitation ratio data. The the-
oretical PDF and CDF are given by (9.5) and (9.7), respectively, with n = 4. Figure
9.2 presents graphical goodness-of-fit analysis: a histogram of the data overlayed
with the theoretical PDF, a comparison of theoretical and empirical CDFs, and a
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Quantile-Quantile (QQ) plot. The graphs show that the fit of the theoretical model
to the data is quite reasonable.

We also examined the assumptions of the model by testing whether the obser-
vations (weekly total precipitation amounts) are distributed exponentially, using the
likelihood ratio test discussed in [14, 19]. The null hypothesis is that the data comes
from an exponential distribution, versus the alternative that the data comes from a
Pareto distribution. Results from the likelihood ratio test indicated that the weekly
precipitation amounts can be assumed to follow an exponential distribution (p-value
= 0.50), with an estimated parameter of ,3 = 0.00648. Figure 9.3 provides his-
togram of the weekly precipitation amounts overlayed with the exponential density
curve with 8 = 0.00648. The graphical fit confirms the results of the goodness-of-fit
test as the density curve closely follows the histogram.
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Additionally, we compared the empirical and theoretical correlation coefficients
between the monthly maximum and the sum. The empirical correlation coefficient
between the sum and maximum precipitation was found to be 7 = 0.871. According
to the results of [20], the theoretical correlation between them should be r = 0.873,
which is rather close to the empirical one.

Our example illustrates that the models involving the ratio of the maximum and
the sum of exponential variables are not only an important theoretically, but are also
promising as stochastic models in actual applications.

Appendix

Proof of Lemma 9. 1. Write the left-hand-side of (9.22) as

(1—g)p ! & j—1 -
T 2 | fenaseeme 02
j=k+2

Note that this is the same as the probability P(T" > k + 2), where T is a negative
binomial random variable given by the PDF

. j—1 a .
s (s+1—1)qs“(1—q)’ GO j=s+ls+2,.

Since P(T > k +2) =1—-P(T <k + 1), we have

00 ] 1 k+1 n 1
s+1 Jj—(s+1) s+1 n—(s+1)
E 1— =1- E 1- )
(s 1 1>q (1-q) (s 1 1>q (1-q)

j=k+2 n=s+1
and the result follows by substituting the right-hand-side above into (9.27). O
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Chapter 10
Least Square Estimation for Regression
Parameters Under Lost Association

Vasudevan Mangalam

Abstract A method is developed to deal with estimating the regression coeffi-
cients when the association among the paired data is partially or completely lost.
Asymptotic properties of the estimators are discussed.

10.1 Introduction

Consider a situation where we have two data sets that are believed to be linearly
related. The independent variable is denoted by x and the dependent variable is
denoted by y. The classical regression analysis gives us least square estimators
for the slope and intercept parameters that are consistent and asymptotically nor-
mal. In this paper the author looks at a situation where the complete data sets for
the independent and dependent variables are available, but the pairwise association
between them is lost. In other words, we have a set of x observations and a set of y
observations without knowing which value of the dependent variable was originally
associated with which value of the independent variable. Such a situation can arise
out of accidental sorting of values that destroyed the association or when the two
data were collected separately or by different people.

The model we work with is the standard linear regression model. Let X,
Xs,..., X, beiid. with mean py and standard deviation ox. Let €1, €2, ..., €,
be i.i.d. with mean zero and standard deviation o, so that €;’s are independent of
X;’s. Finally, let Y; = o + BX; + €;, where « and 8 are unknown parameters. The
observed data consists of {x1, x2,...,x,} and {y1, ¥2, ..., yn} without any infor-
mation on the pairing. In absence of association, it seems natural that we should
define the least square estimator by choosing the permutation of y;’s that would
minimize the error sum of squares. If @(a, ) = mingep, Y 1o (Vri)—(@+pxi))?
where P, represent the set of all permutations of {1,2,...,n}, then our estimators
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of o and § are & and ,3 that satisfy the equation

&, ) = miélcb(a,ﬁ).

10.2 Main Results

In what follows, for any variable x, x,.; represents the ith order statistics of
{X1,x2,...,Xn}

Lemma 10.1. .
(D(&’ ﬂ) = ;IGHPIL Syy(l - rxz;,,(y)) (10.1)

where Syy = Z?:l (yi — )7)2, Ixy indicates the correlation between X and 'y,
and 7 (y) represents the vector obtained by rearranging the values of the vector'y
according to the permutation .

Proof. Let A represent the RHS of (10.1) and let the minimum in (10.1) be attained
by the permutation mp. By the results of standard regression analysis, (see, for
instance, [2]) we know that for any pair of vectors x and y, miny g N [ yi — (a+

ﬂxi)]z = Syy(l—rfy). So for any permutation 7, ming,g Y 1— [V=() — (a—}—,Bx,-)]z
= Sawrm (1 — rf:'r(y)) = Sy(1 - rfn(y))'

4= Sl =riy )

n
= min > oy — (@ + Bxi))?
=1

n
> min min Z(y,,(,-) — (o + Bxi))?
i=1

o,B TeEPy,
= min @(«, B)
a,
= o(@a, p).
Forany 7 € P,,
A < Syy(1— rfﬂ(y))

= min Y iy — (o + Bxi)).

i=1

Thus A < Y7 (Vi) — (@ + Bx;))? Ya and B and Yt € P,. Therefore Ve, f,
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A

A < min > (vre) — (@ + Bx))?
i=1

neP,

= &(a, B).
So A4 < miny g ®(a, B) = P(&, f). O

One of the consequences of Lemma 10.1 is that the permutation that minimizes
@ is not dependent on the values of o and S. If we find the permutation 7z that max-

imizes rf 7(3)” then it minimizes Syy(1 — rf ﬂ(y)) and consequently, ,é = Sxr(y)/ Sxx

and@ = y — BX.

The difficulty is, of course, the computational complexity. If we were to try
out all n! permutations the computational time required explodes at a faster-than-
exponential rate. This problem is tackled by Theorem 10.1. Before that, we need a
preliminary lemma.

Lemma 10.2. Let xy, X3, ..., Xp and y1, V2, ..., Yn be two sets of real numbers
such that x1 < x3 < ..., < Xp, Y1 < V2 < ...,=< Yu. Let P, represent the set of
all permutations of 1,2, ...,n. Then

(1) maxgep, Yty XiVal) = D ey XiVi

(2 minﬂGPn Z:‘l:l XiVr(@) = Z:'lzl XiYn—i+1-

Proof. (1) The proof is by induction. We first prove the result for n = 2.

0 < (x2—x1)(y2—y1) = x1y1 + x2y2 — (X1y2 + x2)1)
= X1y2 + X2y1 = X1)1 + X2)2.

Now assume the result to be true for integers up to and including n — 1. Letting
7 to be an arbitrary element in P,, we will prove that

n n
inyn(i) = inyi-

i=1 i=1
Let j = n(n) and k = 7~ (n) so that w(k) = n. Then

n—1

n
sz'yn(i) = inyn(i) + Xk yn + XnYy;
i=1 i=1
i#k
n—1

= inyn(i) T XkYj ¢t XkYn t Xnyj — Xk Y-
7k
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The expression in the braces, say B, is of the form Z:’:ll X;i yx(;) and hence by
induction hypothesis, B < Zl_l X; yi. Also, applying the result for n = 2 with xy,
Xn, yjand y,, we get X, y; + Xk yn < Xk y; + Xnyn. Hence

n—1

szyn(l) = thyt + Xk Yn + Xny; — XkYj
i=1 i=1
n—1

=Y XiYi + Xy
i=1

n
= Z XiYi.
i=1
(2) Apply Part (1) with y;’s replaced by their negatives. O

Define two perr_nutations Rx and Ry as follows. Ry (i)_is the rank of x; among
{x1,x2,...} and Rx (i) = n — Rx(i) + 1. Let Ry and Ry be defined similarly.
Now define permutations 7r; and m, by m; = R;,IRX and mp = R;,IRX.

Theorem 10.1. Ifif is such that rfn(y) = max{rfm(y), rfnz(y) b then B = Syry)/
Syx and & = y — BX.
Proof. We shall show that

@@, B) = Syy(1 — max{rfm(y), rfnz(y)}),

from which the theorem follows.
From Lemma 10.1, (&, f)=mingep, Syy(1— rfﬂ(y))—Syy(l maxnepn fﬂ(y))

SO it is sufﬁcwnt to prove that maxzep, 7 f a5 = max{ ) s nz (y)} Moreover,

Xﬂl(y
maXgzep, I xyr(y) = max{(maXnEPn rxyr(y)) (mln:rrePn rxr:(y)) }

Note that Z XiVr, () = Z Xn:i Yn:i - Now from Lemma 10.2,
i=1 i=1

neP, n

1 o o
yrrréag; Fxa(y) = |:max - ;(x,-yn(i) —xy):| /sxsy

1 (< _
= - (an:iYn:i —xy> /sty
n i=1
1 n
= (inym(i) —iy'> /SxSy
i=1
= Ixmi(y)-

Similarly, mingep, rxz(y) = Txry(y)- Thus maxzep, r}fn(y) Inax{rm1 )’ mz(y)}.
O
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What we infer from the preceding results is that the least-square estimators are
obtained by ordering the x-values in the ascending order, ordering y-values in
ascending order or descending order depending on which gives a higher coefficient
of determination and finding the standard estimators of the parameters based on this
arrangement. But note that the sign of the slope parameter can be wrongly chosen.
It is possible that a strongly positively correlated bivariate data set yields a higher
coefficient of determination when the variables are ordered in reverse and hence the
method can lead to wrong conclusions on whether § is positive or negative. Thus it
would be prudent to use prior information on the sign of j.

10.3 Almost Sure Limits

In order to find the limits of least square estimators of the regression parameters, we
need the following theorem from [1]. The details of the assumptions are described
in the Appendix.

Theorem 10.2. Let {(Xi(l), Xi(z), e Xi(d)), i=1,2,.. } be a sequence of ran-

domvectors such that foreach j (1 < j <d), {Xl(j), Xz(j), e }forms a sequence

of i.i.d. random variables with continuous distribution function F;. Let ¢ be a real-
valued measurable function on R? such that ¢ satisfies some regularity conditions.
Then

%qu (X,E},?, e X,E‘j’,.)) 2y = E¢ (FT'(U), F5 ' (U), ..., F1(U))

i=1
as n — oo, where U is uniformly distributed over (0, 1).

Note that in the above theorem, no assumption is made about the joint distribution
of X; and Y;. We are now ready to prove the following lemma.

Lemma 10.3. Let F be a continuous distribution function with mean | and stan-
dard deviation 0. Let X1, X2, ..., X, be i.i.d. with distribution function F, and
Y1, Yo, ..., Yy, bei.i.d. with distribution function F. Then,

1 n
_ E an Ynl _)ll.S. //LZ +0,2
n

i=1

and
1
s ,,2 2
; E Xn:i Ynn—it1 —a3 Mmoo —0".
i=1

Proof. Apply Theorem 10.2 with n = 2 and ¢(x, y) = xy, which satisfies the
regularity conditions. Here the limit y is given by
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y=E¢(F'(U). FT'(U))
1
:/ (F~Y(u))*du
0

= /_00 x2dF(x)

o0

=/,L2+02.

The second part of the lemma follows by applying Theorem 10.2 to X and —Y.
O

Lemma 10.4. Let F and G be continuous distribution functions such that there
exists a, b such that G(x) = F(*3%). Assume F has mean p, and standard devi-
ation a1, and G has mean [, and standard deviation 0,. Let X1, X2, ..., X, be
i.i.d. with distribution function F and Y1, Y2, ..., Yy, be ii.d. with distribution
function G. Then,

1 n
n Z Xnii Ynii =4 papa + 0102

i=1

1 n

n Z Xn:i Ynn—iv1 =% pipla — 0107.

i=1
Proof. The result follows from Lemma 10.3 by a linear transformation. O
Next, we look at the asymptotic properties of the estimator. We would hope

that the estimator is consistent and asymptotically normal. Unfortunately, when the
association between x and y is completely lost, we do not get consistency or unbi-

asedness, nor are we able to recover the sign of 8. The limiting behaviour of the
least square estimators are given in the following theorem.

3 a.s. — 2 0_62
Bn =>4 Br =%+ —.
Ox

p =" o =a+ pux (B—P1).

Theorem 10.3.

Proof. From Lemma 10.4,

1
Sxm(y) = ; an:iyn:i — Xy
i=1
—45 E(X)E(Y)+ SD(X)SD(Y)— E(X)E(Y)
= SD(X)SD(Y).

Similarly syz,() =% —SD(X)SD(Y). As 52, =9 V(X) and ,3,, = Sxn(y)/52n
where  is 71 or 7o,
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2 a.s. SD(Y) _ 2 0_62
Pn — so) ~ TVt o
&n = ,)711 - B\nxn
_sa.s. [y — B jix
— at (BB

10.4 Asymptotic Normality

For the asymptotic normality to hold for the least square estimators, we need to make
certain assumptions about the distributions of X and €. In order to make matters
simple, we will not deal here with the case where X and € have general continuous
distributions. We shall assume that both are normally distributed, as a consequence
of which (X, Y') has bivariate normal distribution.

We need to use another theorem from [ 1] as stated below. Once again, details are
given in the Appendix.
Theorem 10.4. Let X; = (Xl.(l), e Xl-(d)) be i.i.d. random vectors. Let F; (1 <
Jj < d) denote the marginal distribution of X l-(j ) \which is assumed to be continuous,
and Fj; (1 < j,k < d) denote the marginal distribution of (Xl-(j), Xl-(k)). If ¢
satisfies some regularity conditions, then

1 n
N p (X;S:Ii)7 S X,Sfll-)) —/n y =4 N0.0% (10.2)
i=1

as n — oo where y is as in Theorem 10.2 and
d 1,y
o? =2 [ [ =@ )dxdy
—170 Jo

1 1
+2 Z /0/0[Gj,k(x,y)—Xy]l/fj(X)l//k(y)dxdy

1<j<k<d

where G jx(x,y) = Fjx (Fj_l(x), Fk_l(y)).
Lemma 10.5. Let (X1,Y1), (X2,Y2), ..., (Xu, Yy) be i.i.d. bivariate normal with
each component being N (0, 1). Then,
1. \/L; Z?:l (Xn:i — Yn:i)2 =70
2. =Y [XniYni = Xn¥u — 1] >4 N (0,72%)
where 12 = % [E (XZYZ) + 1].
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Proof. For Part 1, apply Theorem 10.4 with n = 2, ¢(x, y) = (x — y)2. It can be
verified that when the distributions are normal, the conditions of Theorem 10.2 and
Theorem 10.4 are satisfied. It is easily seen that for this case, the asymptotic mean
and variance are both zero, and hence the result holds. As for Part 2,

2Xn:i Yni = X,%;,' + Ynz;i - (Xn:i - Yn:i)2 s

SO

n n n n

2X:Xn:iyn:i = ZX,Z + Z Y,'Z - Z(Xn:i - n:i)2

i=1 i=1 i=1 i=1
1 < - — 1 &[X2+7Y2 — —
WE[Xn:iYn:i_XnYn_l]z%;[%_1]4_\/5)("}/"4_017(1)

from Part 1. By the Lindeberg-Lévy central limit theorem (See [3], for instance),
the first term of RHS goes to normal with mean zero and variance equaling

X2 +v?
()
2

[V (X?)+V (Y?) 4200V (X?,Y?)]

N — ] — A —

[24 cov (x>, Y?)]

[E(X?Y?) +1]

N

)

ﬁ X, Y, —P 0because /1 X, is asymptotically normal, and hence O (1), and
Y, =2 0. The result now follows from Slutsky’s theorem. O

2
Note: The asymptotic variance 2 is equal to V(f ) if X;’s are independent of Y;’s,
whereas t2 =V (Xz) if X; =Y, foralli.

Lemma 10.6. Let (X1,Y1), (X2,Y2), ..., (X,,Yy) beiid. bivariate normal such
that X1, X2, ..., Xy areiid N(uy,01)andYy, Ya, ..., Yy beiid N(uz,02).
Then,

n
% Z [Xn:i Yoi — (Yn?n + 0102)] _>d N (07 772)

i=1
2.2 2 2
where n? = 01202 E |:(X;1‘“) (Y;é“) + 1] )

Proof. LetZ; = Xizl and W, = Y’U;z“z By Lemma 10.5,

a1

1 <& o
%Z [Zn:i Wn:i — Zan — 1] —)d N (O, n2t) )
i=1
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The left hand side is equal to

n J—
«/ﬁ;wz i; [X,,;,-Yn;,- - (YnYn + 0102)] and hence

12022 [E (ZZ W2)
+1] which equals 7. |

We are now ready to prove the asymptotic normalities of ﬁn and @&,.

Theorem 10.5. If (X1,Y1), (X2,Y2), ..., (X,,Yy) are ii.d. bivariate normal,
then the least square estimators of the regression parameters under lost association
are asymptotically normal.

1. Vi (B = 1) =7 NO.)
2. Jn(dy —ay) =4 N(O,")

where By and oy are as in Theorem 10.3

Proof. Bn = sx,,(y)/s)%n where 7 is m; or . We shall prove the case of 7 = 7.
The other case is similar. (It may fluctuate between the two permutations giving
positive or negative values for ;. In that case the subsequences will converge to 81

or —f1). As 52, =45 o2,

i (B = 1) = i (B2 - 202

2 2
Sxn Ox

1 ¢ -
=n (; > XwiYmii — xy/s;‘in)

i=1

J_UZZ Xn:i Yo — (_Yn+0102)]
X

i=1

The asymptotic normality of ,3,, now follows from Lemma 10.6. The asymptotic
normality of &, follows from that of ﬂn and the factthat & = y — ﬁx |

10.5 Partial Loss of Association

It is conceivable that only for part of the data the association between x and y is
lost. For the rest, we may have the full data set with the one to one correspondence
intact. In such situations we expect consistency and asymptotic normality for the
least square estimator to hold provided the proportion of observations for which
association is lost tends to zero.

As this is a rather severe restriction, the author suggests alternative estimators
for B and « as follows. Let s2, be the standard estimator of the error variance, s2,,
be the standard estimator of the variance of the x variable, and 8 be the standard
estimator of 8. These estimators are calculated using the part of the data for which
association is not lost.
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- R 2
B = sen(y)[Bn” — .

Assuming that the number of observations for which association is intact tends
to infinity as the sample size tends to infinity, this new estimator is consistent. This
is a straight forward consequence of the previous results. Its asymptotic normality
is, at present, not as clear.

10.6 Conclusion

There is scope for further studies on this topic. Investigating MLE’s is one possible
extension. Another is to generalize the idea of lost association. In general, loss of
association does not have to be a total hit or total miss. It is possible that for each
Xx;, we have a set of y;’s that x; may be associated with, and this set may range
from a singleton to the entire set of y;’s. On one extreme we have full association
information and on the other we have none. Least square estimation and maximum
likelihood estimation for such a general case can be a subject of future investigation.

Appendix

The content of this appendix is an excerpt from [1].

Let {(Xi(l), Xi(z), e, Xl-(d)), i =1,2,...} be asequence of random vectors such
that for each j (1 < j < d), {Xl(j), Xz(j), ..., forms a sequence of independent
and identically distributed (i.i.d.) random variables with distribution function Fj.
Let X,Ej-) denote the i th order statistic of {Xl(j), Xz(j), ... X,Ej)}. Define, for0 < ¢ <

N

1, F~(¢t) = inf{x : F(x) > t} for a distribution function F.

Let ¢ be a real-valued measurable function on R Letx = (x1,...,xq) for
0<x,x1,...,xq < 1.Define
Y(x) =¢ (F{ '(x1)..... F; ' (xa)), (10.3)
y(x) = ¥(x,x,...,x), (10.4)
oY (x i

vi(x) = #Rx ,,,,, X, 1=<j=<d (10.5)
Xj
0%y (x )

Vjk(X) = 3 id ), <jk<d. (10.6)
X j 0Xk

We introduce conditions on ¢.
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(C1) The function v (u) is continuous atu = (u,...,u),0 < u < 1. Thatis, ¥ is
continuous at each point on the diagonal of (0, 1)¢.
(C2) There exist K and c¢ > 0 such that for (x1,...,x4) € (0, co)d U (1—=co, l)d,

d
W, xa)l < K[ 1+ Iv(x))]

Jj=1

(C3) The functions, [x(1 — x)]3/2[1p,- X2 (1 < j < d),and [x(1 — x)]?/?
[¥jk(x,...,x)| (1 < j,k <d)are Riemann integrable over (0, 1).
(C4) For all large m, there exist K = K(m) > 1 and § > 0 such that

[ (y) =¥ () =y —x. VY (x))|
d
<K Y10 =00k =) 1+ ¥l

J.k=1

ifx = (x,....x),y = (V1,....y4) € (0,14, [ly — x||¢, < &, and for
l<j=d yj(d=y;)>x(1 —x)/m. Here|yl¢ := |y1] + -+ [yal
denotes the £;-norm of y and V/(x) the gradient of .

Theorem 10.6. Let {(Xl-(l), Xl-(z), e, Xl-(d)), i = 1,2,...} be a sequence of ran-

dom vectors such that foreach j (1 < j <d), {Xl(j), Xz(j), ..., } forms a sequence
of i.i.d. random variables with continuous distribution function F;. Suppose that
¢ satisfies the conditions (C1)—(C2), and that y, as defined in (10.4), is Riemann
integrable, then

1 " ) @)\ a-s. -
;Z(p(Xn:i""’Xn:i)—_) 7
i=1
asn — oco. Here j = [y y(y)dy = E¢ (F{'(U), F; ' (U), ..., F; (U)) and
U is uniformly distributed over (0, 1).

Theorem 10.7. Let X; = (Xl.(l), e Xl-(d)) be i.i.d. random vectors. Let F; (1 <
Jj < d) denote the marginal distribution of X l-(J ) which is assumed to be continuous,

and Fjr, (1 < j, k < d) the marginal distribution of (Xl-(j), Xl-(k)). If ¢ satisfies
conditions (C1)-(C4), and that y, as defined in (10.4), is Riemann integrable, then

1 é _ dis
NG Y ¢ (X,E;l,-)’ s Xﬁfl,-)) —Vn7 5 N, 6% (10.7)

i=1

as n — oo where y is defined as in Theorem 10.6 and
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d 1 y
02 =23 /0 /0 * (1= ) ()W ()dxdy
j=1

1ol
+2 Z /0/0[Gj,k(X,Y)—Xy]l//j(x)l/fk(y)dxdy

1<j<k<d

where Gj(x,y) = Fjx (Fj_l(x), Fk_l(y)).
If we arrange the random variables in a tabular form as below:

Xl(l) Xl(j) de)
Xz(l) sz Xéd)

In Theorem 10.6, we just require i.i.d. for each column and no further assumptions
are made on how the columns are related. We need a stronger assumption in Theo-
rem 10.7, namely, the rows are i.i.d. random vectors. Interestingly, the variance of
the limiting normal only depends on the 2-dimensional marginal distributions.

We collect some remarks about the various conditions on the function ¢.

Remark. (i) Conditions (C1) and (C2), in general, are easy to verify.
A sufficient condition for condition (C4) to hold is that the second par-
tial derivatives of ¥ are continuous, and there exist K and @« > 0 such

that [Y 4 (y1.....ya)| < K X0y Wk (... yo)l and [k (x. ... x)| =
O(x(1—=x)]7%) for1 < j,k <dasx — 0+ orasx —> 1—.

(i) By a compactness argument, Condition (C1) is shown to be equivalent to

(C1”) For any ¢ € (0, %) limg_,q w(c,8) = 0 where

w(c,8) == sup|¥(x1,...,xq) —y(¥)| (10.8)

and the supremum is taken over all ¢ < x;,x2,...x4,y < 1 — ¢ such that
|xi —y| <38.

(iii) Let ¥(x,y) = [(x +y)/2]*[1 — (x + y)/2]%. 1t is easy to verify that for
a > —1/4,  satisfies conditions (C3).
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Chapter 11
On Tests of Fit Based on Grouped Data

Sherzod M. Mirakhmedov and Saidbek S. Mirakhmedov

Abstract The problem of testing the goodness-of-fit of a continuous distribution
for a set of n observations grouped into N equal probability intervals is considered.
It is assumed that N — oo as n — oo. Let 1, ...,ny be the numbers of obser-
vations in the intervals. We show that within the class of tests based on statistics of
the form f (n1) + --- 4+ f (nn) the classical chi-square test is optimal in terms of
the Pitman’s and the Kalenberg’s intermediate asymptotic efficiencies but it is much
inferior to tests satisfying Cramer condition in terms of the Kalenberg’s strong inter-
mediate and the Bahadur’s exact asymptotic efficiencies. For the chi-square statistic,
a probability of large deviation result, likely to be of its own interest, is proved.

11.1 Introduction

The classical goodness-of-fit problem of statistical inference is to test whether a
sample has come from a given population. Specifically, we consider the problem
of testing the goodness of fit of a continuous distribution F to a set of n observa-
tions grouped into N equal probability intervals. A large class of the tests based on
statistics are of the general form

N
Ry =" f(no. (11.1)
k=1
where 11, ..., ny are the numbers of observations in the intervals. In the sequel the

statistics R{, is called f-statistics, a test based on f-statistics is called f-tests. Three
most popular cases of f-statistics are
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N
2 2
Xy =2
k=1

is called the chi-square statistic,

N

Ay =) mlnm
k=1

is called the likelihood ratio statistic,

N
tr =Y A inm=r}
m=1

is a number of intervals containing exactly r observations, here f} {A} is the
indicator of the event A. Particularly p¢ is known empty boxes statistic (see, for
instance, [16]).

We consider the problem of testing the null hypothesis Hy: f(x) = F'(x) = 1,
0 < x < 1, versus the sequence of alternatives

Hy: f(x) =1 +d8n)l(x), (11.2)

where constant d > 0, [(x) is a known continuous function on [0,1] such that

1 1 1
/ I(x)dx = 0, / I?(x)dx =1, and / [1(x)|? dx < oo,
0 0 0

8(n) — § < oo will be chosen so that the power for a f-test of size w has a limit in
(w,1). The problem of testing Hy against H; is called problem (Hy, Hy).

Holst [4], Ivchenko and Medvedev [6], and Gvanceladze and Chibisov [3] have
shown that for the problem (Ho, H,), for §(n) = n~/2 the power of the f-tests
tends to the significance level as n — oo whenever N — oo; hence such alterna-
tives can not be detected by f-tests. In [20], see also [30], it was pointed out that if
we let N — oo then a sequence of alternatives that convergence to uniform must
be in the form (11.2) with 8(n) = (na)~'/*, where @ = n/N, in order to keep
the power bounded away from the significance level and unity, hence the f-tests
do not discriminate alternatives (11.2) with §(n) = o ((n)~'/#). This is poor in
comparison with other tests based on empirical distribution functions, for example
the Kolmogorov—Smirnov and Cramer—von Mises tests, which can detect similar
alternatives at distance O (n_l/ 2). On the other hand not always we need to con-
sider the alternatives converging to hypothesis with the extreme rate of O (n_l/ 2).
Moreover, concerning the choice of the number of groups in chi-square test there is
a well-known result by [17] stating that the optimal number N increase with n as
N=0 (nz/ 5 ) Hence it is unnatural to keep fixed number of groups when number
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of observations goes to infinity. Nevertheless, because of §(n) = (na)~'/* the
number of groups should go to infinity slowly in order to discriminate alternatives
converging to the hypothesis with the rate close to O (n~'/2).

We are concerned with asymptotic results when N = N(n) — oo as n — oo.
This case has been studied intensively by many authors. We refer to [1, 4, 6, 9—

11,15, 18-20, 27, 29, 30]. For Rf, [7,8,21,36] prove: the central limit theorem
under mild condition together with Berry—Esseen bound, Edgeworth type asymp-
totic expansion with exact formula for the first three terms, specified for statistics
X 12\,, Apn and p,, and Cramer type large deviation result under Cramer condition
Eexp{H |f(§)|} < oo, IH > 0, where £ is the Poisson(«) r.v. with @ = n/N.
We refer also to [34] where under Cramer condition the Chernoff type large devi-

ation result for R 1{, was proved. Note that the statistics Ay and p, satisfy Cramer
condition whereas the chi-square statistic X 1%, does not. Nevertheless [30] obtain
asymptotic results for Chernoff type large deviation probabilities of X 1%, and Ay.
These probabilistic results have been used to study asymptotic efficiencies (AE) of
the f-tests. Details of the corresponding results are as follows.

There are two basic ways of comparison of tests. One of them is in principle
based on asymptotic analysis of the power of the tests. A test having maximal
power within a class of tests under consideration is called asymptotic most pow-
erful (AMP) test. AMP test is also first order efficient test, and it may be not unique.
In such a case an asymptotic behavior, as n — 00, of the difference in powers of two
AMP tests is of interest; this situation gives rise to the concepts of second order effi-
cient tests. The definition of the second order efficient test adapted to our problem
is given later.

Another method of comparison of two tests of the same level is based on com-
parison of the number of observations needed to get same asymptotic power, when
number of observations increases. If we have two tests with corresponding numbers
of observations n; and n,, then the limit of ratio n,/n; is called the asymptotic
relative efficiency (ARE) of test 1 with respect to (w.r.t.) test 2. To investigate AE
of a test we consider that ratio where n; and n, corresponds to that test and the
AMP test respectively. AE of a test depends on three parameters: the level w,, the
power f3,, and the alternative H;, which may depend on n. When letting n go to infin-
ity, three concepts arise: Pitman approach when w, — @ > 0, H; — Hj in such
rate that 8, — B € (w, 1); Bahadur approach when v, — 0, 8, — B € (0,1), H;
is fixed, i.e., does not approach the hypothesis; Kallenberg intermediate approach
when w, — 0, 8, — B € (0,1), Hy — Hy but more slowly than in Pitman
case. Optimality of a test can be expressed by first order efficiency, which means
that ny(w, B, H1)/n1(w, B, Hy) converges to 1, where the limit is taken accord-
ing to the efficiency concept involved. The problem of finding of the limit of ratio
na(w, B, Hy)/ni(w, B, Hy) is a very difficult problem as it usually reduces to find-
ing of the ratio of what is called slopes of the tests under consideration, see, for
instance, [5, 28].

Let £ be the Poisson(a) r.v. with @« = n/N; P;, E;, Var; stands for the proba-
bility, expectation and variance under H;; A; y and ol% ystands for the asymptotic
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value of E,-RI{I and Var,-Rj{,, i =0, 1, respectively. Put
gE) = fE —EfE —yE—a).y =a cov(f(§).§) (11.3)
o*(f) = Varg(§) = Var f(§) (1 —cor® (f(§).§)) .

From Theorem 2 of [21] it follows that if

_ Elg®

= 28N o, 11.4
KN NG - (11.4)

asn and N — oo, then

7 1
Vi = sup| Py {Rf < xouy + Aiw | — 9()] = O (KN + W)’ (11.5)

where @(x) is the standard normal distribution function.

Remark 11.1. The statistics Xj%, and Ay satisfy the condition (11.4) if and only
if na — oo. But for the statistic u, (11.4) is valid under additional conditions
for r and o, namely, if « — 0 and no — oo then (11.4) satisfies for u, with
0 < r < 2;if « is far away from zero then (11.4) is still true for u,, r > 0, if
o« —InN —rInlnN — oo.

In what follows we will assume that noe — oo.
Asymptotically Most Powerful Test. It is known, see [4,7,8], that under alternative
(11.2) with §(n) — 0

Aoy = NEf(£),02y = No2(f),0ly = oay(1 +0(1)), (11.6)
v ()L Ain(f) = Aow (f)) /VNo(f) = \/?SZ(n)p(.ﬁ a)d? (1+o0(1)),

(11.7)
with p(f &) = corr (f(§) — y§.6% — 2o + D).

Let S(f) be an asymptotic power of the f-test of a size w. If (11.4) is satisfied
then (11.5) and (11.7) imply

B(f) =@ (@Sz(n)p(ﬁ a)d? — uw) iy = P71 —w). (11.8)

Hence the functional p( f, @) plays a key rule in determining of the asymptotic
quality of the f-test. Its meaning is clarified by the following (see Lemma 1 by [8])

o(f. @) = lim corro (R,{,,X},). (11.9)
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Hence |p(f,@)] < 1, and |p(f, )] = 1 for any « only for chi-square test. The
equality (11.8) means that f-test does not detect alternatives (11.2) with §(n) =
o ((na)=14).

In (11.2), let §(n) = (na)~/*. Then for the problem (Ho, H,) the chi-square
test is AMP within class of f-tests for any «. Nevertheless if @ — 0 or « — oo then
there exist other AMP tests also, because in these cases we may have p( f, o) — 1.
For example, if o« — 0 and A? £(0) # 0, where operator Af(x) = f(x+1)— f(x),

then s
~ 22O 6 02);

p(fia) =1 6 A27(0)

if & — oo for the statistic Ay

p(fie) = 1= o (1+0(1).
o

Second Order Asymptotic Efficiency. Comparison of the AMP tests, when o — 0
or ¢ — 00, can be based on the notion of the second order efficiency. Set =
d?/v2 — ug,, where w is a size of f-test and ug, from (11.8), By (R{,; 1) stands
for the power of the f-test of a size w. Ivchenko and Mirakhmedov [7] show the
following asymptotic expansion of the power 8, (X 1%,; ) of chi-square test

Bu(X3:0) = @ () + en(X3:9) (1 4+ 0(1)),

where

exp {—92/2} (1 - 92 9d? ne n
W(X3:0) = +—+ V25 (? —+—)),
i) = e 3y T2 UV T2

ifa — 0,

2 2
ey = SO (0
27N V2
if ¢ — oo.

Here S;(x) = —{x} 4+ 0.5, {x} denotes the fractional part of x. The func-
tion S1(x) is well known in the theory of asymptotic expansion of the cumulative
distribution function of lattice random variables (r.v.’s). It arises here because the
chi-square statistic X 1%, is a lattice random variable with span equal to 2.

Definition. The AMP f-test is called second order asymptotic efficient (SOAE)
with respect to chi-square test, if its power has the asymptotic expansion

Bu(RY.9) = @ (9) + ea(f39) (1 + 0(1)),

with g,(f: ) — 0and &,(f: ) = ex(X3%: ) (1 + o(1)).
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The conclusion of [7] is as follows. Let « — 0, then there exist SOAE f-tests
only if « = O ((na)~Y/2),i.e., n = O (N3/*). For example empty cells test based
on (o is SOAE, but likelihood ratio test is not SOAE. If « — oo, then SOAE does
not exist.

Pitman Efficiency. Under certain regularity conditions (see, for example, [2]), the
efficacy of a test based on statistic, say V, is given by e(V) = u%, / 0%,. Here py and
0%, are the mean and variance of the limiting normal distribution under the sequence
of alternatives when the test statistics has been normalized to have limiting standard
normal distribution under the hypothesis. In such a situation, the Pitman’s ARE of
one test with respect to another is the ratio of their efficacies.

Because of (11.6) and (11.8) the alternatives (11.2) with §(n) = (ne)~"/* are
form P,;-the family of Pitman alternatives: f-test of a size w,(f) — @ > 0 and
p(f, a) is far away from zero has the power (see (1.8))

Bu(f) = B() = & (272d%0(fe) — o) € (@, 1).
For the efficacy e(f) of the f-test we have

e(f) = x5 (f) =271d**(f ).

Hence the Pitman asymptotic efficiency of f-test is determined by functional
p(f, ) and within class of f-tests the chi-square test is asymptotic most efficient
(AME) in Pitman sense; the Pitman efficiency of f-tests goes down as number of
intervals N increases for a given sample size n. These results have been proved by
[4,6,20,30].

Bahadur Efficiency. Another extreme family of alternatives is B,,-Bahadur (as
well as Hodges—Lehman) family of alternatives when alternatives do not approach
the hypothesis, i.e., §(n) is constant. The Bahadur’s AE of f-tests in the family
B,i; have been developed by Ronzhin (1984) who showed, for a certain subclass
of f-tests, that whenever B, (f) — B(f) < 1, —n~'log w,(f) converges to limit
(which is called slope of the f-test to the alternative) of specifies the Bahadur AE of
f-test. This limit is determined by the logarithmic rate deviations probabilities (for
deviation of order O(+/N)) under Hy, which require restrictive Cramer’s condi-
tion (see (11) below) on the test statistics. In particular, this condition excludes the
chi-square statistic. A comparative analysis of chi-square test’s Bahadur efficiency
relative to the likelihood ratio test was carried out by [30]. They showed that likeli-
hood ratio test is much more Bahadur efficient than the chi-square test, in contrast
to their relative Pitman efficiency. In a similar setup, the ARE, as « — oo, of the
chi-square and likelihood ratio tests were studied by [15] based on the analysis of
probabilities of large deviations of various orders.

Kallenberg Intermediate Efficiency. The situation when §(n) — 0 but slower
than that in the P, give rise to the Kallenberg’s intermediate family of alternatives
of three types: K-family of alternatives (11.2) with §(n) — 0, §(n)(na)'/* — oo;
K ¢-subfamily of K,; with 8§(n) = o ((naz)_l/G) if o > 1, and §(n) =
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0 ((na)_1/6) ifa < 1; K g -subfamily of K,y with §(n)=0 (not)_l/4 logl/4 N).
Actually such division of family of intermediate alternatives becomes from prob-
ability of large deviations results presented below in Sect. 11.3 and because of
relation (11.7). Following the logic of Bahadur’s approach, intermediate AE (IAE)
(between Pitman and Bahadur settings) of f-tests for the family of intermediate alter-
natives (Ku, K176 and K J@) can be measured by the logarithmic rate of decrease
of the test size when the power is fixed. Therefore, [8] consider as a measure of the
performance of f-test, the asymptotic value of a slope

¢®(RL) = —log P, {R{, zNAlN(f)}. (11.10)

ARE of one test relative to another is defined as the ratio of its asymptotic slopes.
For Pitman’s alternatives this is equal to the Pitman’s asymptotic relative efficiency,
whereas for intermediate alternatives it is related to the IARE in weak sense intro-
duced by [5]. Ivchenko and Mirakhmedov [8] extended this above said result on the
Pitman’s and the Bahadur’s efficiencies properties of the chi-square test: chi-square
test is still optimal within the class of f-tests in the Kallenberg’s weak IAE (i.e., for
the family K /5;) but in the Kallenberg’s strong intermediate efficiency (i.e., in the
family K,y except Kj/e) it is much inferior to those statistics satisfying the Cramer
condition, particularly to likelihood ratio test.

It follows from what has been said above, that the chi-square test is AMP, SOAE,
and AME in the Pitman’s and the Kallenberg’s weak IAE senses, but it losses opti-
mality property in terms of the Bahadur’s and the Kallenberg’s strong efficiency. For
the f-tests satisfying Cramer condition AE for all range of alternatives (11.2), i.e.,
for family of alternatives P,;, K,; and B, in the situation when « is far away from
zero and infinity have been studied also. AE of the chi-square test in the family of
alternatives K/ has been an open problem.

Our main purpose here is 2-fold. On the one hand we wish to cover that existing
gap in the study of AE of the chi-square test; with this aim we prove a probability
of large deviation result, likely to be of its own interest. On the other hand we will
extend the result of [8] regarding to AE of chi-square test in the family K,;; except
K¢, for the cases « — 0 and o — oo.

11.2 Main Result

We assume na — o0. Also we continue to use the notations as in (11.3), (11.6),
(11.7) and (11.10). The main conclusion of the present paper is the following.

Theorem 11.1. /. If0 <co <o <c¢1 < o0 and

Eexp{H | f(§)|} < oo, (11.11)

for some H > 0, then in the family of alternatives K .
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w(R ) d4

iy = g P U@+ o(),

2. In the family of alternatives K¢

w(X ) d4
P = (o).

3. In the family of alternatives Ky if

83(n)(na)?log™' N — oo, (11.12)
then o(x2)
ey (X .
nadt(n) o).

This Theorem 11.1 together with the results of [7, 8, 30] implies that chi-square
test is optimal within class of f-tests in Pitman AE and Kallenberg IAE senses but
it is much inferior to those statistics satisfying the Cramer condition (particularly to
likelihood ratio test and w,,r > 0, tests) in Kallenberg strong IAE under condition
(11.12) and Bahadur AE senses. These covers almost all range of alternatives. The
only remaining gap in the study of the intermediate efficiency of the chi-square test
is in the family K,; with

c2(na?)™V8 < §(n) < c3(na)™V0log"? N

fora > 1,
and

cz(not)_l/6 <én) < C3(n0l)_1/6 logl/3 N
fora < 1.

Remark 11.2. An alternative approach to testing of uniformity [0,1] is to construct

tests based on “spacings”. Let X1, < X2, < ... < X, , be the order statistics
of the sample X1, Xa..... Xp, Wb = Xpsn — Xm—tys»m = 1,2,....N’,
W(s) = 1 — Xnvs,4, with notation X, , = 0 and X,41,, = 1, be their s-

spacmgs N =[mn+1)/s], N=N"if (n+1)/sisanintegerand N = N’ + 1
otherwise; W = (Wl(sn), e,
with n, but s = o(n).

WA(,SL) The step of the spacings s may increase together

The order statistics Xon, X1n, ..., Xn,n, Xn+1,n divide interval [0,1] to s + 1
subintervals (groups), that is we again, actually, deal with method of grouping data.
In contrast to above considered method here the ends of intervals are random and we
are using, for a statistical procedure, the length of intervals instead of frequencies of
intervals. Most common among tests based on spacings are tests based on statistics
of the form
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N
Rewy =3 1 (0 +1w8)).
m=1

AE properties here alike to those of f-tests (the step of spacings s plays the role
of «). For example: such tests can detect alternatives (11.2) with §(n) = (ns)~¢,
e € (0,1/4], AMP test for the alternatives (11.2) with §(n) = (ns)~'/4 is the
Greenwood’s test based on statistic

N
G =Y (n+ 1)W,§j3)2.
k=1

While considerable attention has been devoted in literature to type R 1{, (W) statis-
tics, we are not in position to give here all the details of existing results. Reader can
find detailed information, applications, and references, for example, in papers by
[11,13,23-25,32,33,37]. We wish only to refer to [9—11] where the Pitman’s ARE
of chi-square test and Greenwood test with s = [«] were studied. They have shown
that if s — oo then these two tests have same Pitman efficiency, but for fixed s (that
corresponds to the case 1| < a < ¢ < 00) spacings tests are preferable to compara-
ble chi-square procedures. From Theorem 2.1 and results of [26] it follows that the
same results are still true for Kallenberg IAE of chi-square test and spacings based
tests.

Proof of Theorem 11.1 For proof of Part 1, we refer to [8]. To prove of Part 2 we
note that

e?(RY)

—log Py {RI{, > xn(f)o(f)VN + Nf(é)}

f_
—log Py { Ry —NJG©) | g82(n)p(f,a)d2(l +0(1))}

o(/INN

because of (11.10). Therefore, Part 2 follows from Theorem 11.1 of Sect. 11.3,
using the facts that p( f, @) = 1 for the chi-square statistic and that — log @(—x) =
271x2(1 + 0(1)), as x — oo. O

Proof of Part 3. We use an approach similar to that of [30]. We will write n ~
B(k, p) to mean that a r.v. 5 has the Binomial distribution with parameters n and p,
0<p<l;andv(n) = [a + Vo + dznot(gz(n)] + 1 where [a] is an integer part
of a. We have

P{X3 >NA) =P {XJ%, —n@+1)> xN(_f)vzna}

> ((hm — @) — @) > d*nas*(n) (1+0(1))§

N
m=1
N

> (O — @) —a) 20/771 = v(n)

m=2

=P
=P P {m =v(n)}
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N—-1
=P ((im—a)*—a) 20} Pin=v)}.  (11.13)
m=1

Here 7, ~ Bi (n —v(n), (N — 1)_1). Puta = (n —v(n))/(N — 1). It easy to see
thatv(n))/n = (N7' + dS(n)N~1/2) (1 + o(1)) and @ = a(1 + O(N ! +dé(n)
N~1/2)). We have

N-1 N—-1
PIY ((m—)?—a) = 0} > P> (im—&)? = (N - 1)a}
m=1 m=1
N-1
=P ((lm—@)?*—a) = (v(n) —a)
m=1
Yot (Gim —8)2 = &)
=P ds 1
V26— - e
> ¢ >0, (11.14)
because of (n — v(n))a = no (1 +o0(l)) — oo, and hence the Central Limit

Theorem for chi-square statistic ZZ;% (fm — @)? is valid, see above Remark 11.1.

Set g(x,p) = xlog(x/p) + (1 — x)log((1—x)/(1 —p)), x € (0,1) and
p € (0,1). The following Lemma 11.1 is quoted from [30]. O

Lemma 11.1. If§ ~ Bi (k, p) then for integer kx
P& =kx} > 0.8 2rkx(1 —x)) Y2 exp{—kg(x, p)}.

Note that 1 ~ B(n, N~1). Therefore due to Lemma 11.1 we have

P{nm=vin)}=>c [v(n) (1 — v(n)n_l)]_l/2 exp{ —v(n)log(a™'v(n))

—n(1 —n"v(n))log %}

> ¢ (v(n)) "% exp {—v(n) log(a@™ v(n))} .
Hence
_log P im =v(m)i _ clog v(n) +v(n)log(a~"v(n))
nad4(n) - nos*(n)

a+8(n)/no . v(n)
c——————log —

nad*(n)

1 1
=¢ |:7’l84(n) + 83(’1)\/@] log N =o0(1), (11.15)

because of (11.12). The Part 3 follows from (11.13),(11.14) and (11.15).
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11.3 Large Deviation Results

We recall from the notations of Sect. 11.2: £ is Poisson () r.v., « = n/N,y =

o~ leov (f(§).6),

g&) = fE& —EfE) —yE—a) .o (f) = Varg(§)
= Varf(€) (1 —corr* (f(£),£)). (11.16)

The following result on the large deviation probability of the chi-square statistic
stated in Theorem 11.2, is likely to be of its own interest.

Theorem 11.2. Ifa > 1 thenforallx > O0and x = o ((n/ot)l/ﬁ), if o < 1 then for
allx >0andx = o ((not)l/ﬁ) one has

P {XJ%, > xv/2na + n(o + 1)} — (1—®(x) (1 +o(1)).

Proof. Let &1, &, ... be independent r.v.’s with common Poisson («) distribution.
Also let Cg (§) be a cumulant of the kth order of ar.v. £ and

SN—Z(sm @), XIN—Z(nm— X%N—Z@m—a) 0

Lemma 11.2. For any fixed k > 2 and large enough n
Ce(X7 y) = Ce(X7 ) (1 + 0 (1))

Proof. 1t is well known that L((i11,...,0n8)) = L((&1,....EN)/SN = 0),
where £(X) denotes the distribution of the random vector X . Hence

E(Xy)F = E ((X2)"/Sy =0). (11.17)
On the other hand E ((Xlz,N)ke”SN) =E {ei’SNE ((XIZ’N)k/SN)}. Integrating

w.r.t. T both side of this equality over interval [—m, 7], and taking into account
(11.17) we have

k Tk Sy
E (X? =d E (X? exp{ir—} dr,
( l,N) n —_ ( l,N) «/ﬁ
where
1 nle"

dy = (2m/nP{Sy =0}) " = P
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Hence, putting §m = §,, — a we have

E(X2 )k d Zii[”ﬁE[gk‘ §2klex {irSN}]dr

o n1=1 K 1 Jmn At O Vn ’
(11.18)
where Y} is the summation over all I-tuples (k1, ..., k;) with positive integer com-
ponents such that k1 + --- + k; = k; Z;’ is the summation over all
I-tuples (J1, ..., j;) with components not equal of each others and such that j,, =

L2,...,Nym=1,2,...,1
Putting Sy; = Zzl‘=1 &, we have

wn B B S
2k 2k . N
/_nﬁE[Eh‘-...-Sjllexp{ztﬁ”df
T . N Sn—S
_ gleza . g { N,IH[E { N N,l}
/;nﬁ |:E“ é]/ exp lr—ﬁ exp lr—ﬁ
2 -1 w/n 2 -1
_ (1—L) d7:+/ exp - (I—L)
2 N - 2 N

<[ (owfie <) Jare £ 8]

— exp

w/n 2 i -1
x/ expl - o (1 - —) ™ 5 1+ U5, (11.19)
./ 2 N
We have
g23 2
E exp l:/gg’} ‘ =exp{—2asin2 Zj/ﬁ} §exp{—%r2} , (11.20)

since sin? u/2 > u?/n?, |u| < 7. Put A, = 3N~Y2 4 (na)~1/2, then /nVar3/?
Em/E |Em|® = A, !, Taking into account this inequality and using Assertion 11.4
and (11.20) by some algebra we have

g z . SN — SNy
< E[8R. gk {/ E { —}
= [E“ i ] ezeant | L

72 I\t
_exp§—7(1_ﬁ) } ar
Sy —S
+/ (‘Eexp{it—N N’IH
7w /nzlt]2(64,) NG
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? 1\ k g2k
+exp{—7(l—ﬁ) dty < C4AL E [Szllu..-*;‘jll].
(11.21)
Lemma 11.3. For any integer v > 2 one has
B lv/2]
EE, =) cdl (11.22)
with
O0<e,<1,1=12,...,[v/2]. (11.23)

Proof. We have Ee'™ = exp {o (¢'® — 1)}. Applying here the Bruno’s formula
we find

v [v/2]
- 1
Vol ko+...+ky
EE V.ZO( U '(m')km =! ch Voz
m=2
where ) is summation over all non-negative k», . . ., k, such that 2k, +. ..+ vk, =

vand! =k, + ...+ ky,cip =Y [Lnhes W Lemma 11.3 follows.

In particular, from Lemma 11.3 it follows that EEX*+1 < CEE? and EE2k+2 <
Cok2aEE?* . Using this fact after some algebra we obtain

1 2k 22k; @2 0 2 72 ) !
|J2|<—E[§ 1'---'§j1151v,1]/ Thexp) —— 1_N dt

—00
ak?l + k212 [~ .
<C———E [Zj.l"js Zjl"g] (11.24)
It is obvious that
I
-2 E[Zkl-... 52"’](1+0(N)). (11.25)

Apply (11.21), (11.24) and (11.25) in (11.19) to get

[ (e et e

2 3
=@(1+0(k7+%+%+\/#_))12[52’” -...-éj.l"l]. (11.26)

Using Stirling’s formula it is easy to see that d, = (27[)_1/2 (1 + O(n_l)).
Inserting this and (11.26) into relation (11.18) we have
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3
E(X3y) = E(X2y) (1 +0 (% + ﬁ + J%)) .12

Lemma 11.2 follows from Assertion 11.3, see Appendix, and (11.27), because
Ce(X3) = Ce(XT y) -
Let @ > 1. Due to Lemma 11.3 and Stirling’s formula we have: for k > 3

£ (6282

k—2 ~ _ ~
< (k)2 23+ ok < (k1) (21°k1/<k—2>a) Varg2 < (k)2 (2'%a)* > Varf2.

k
= zkEgr%,k < (2k)12* Zal < (kN2 2L 2¢ (Zk)
I=1 (k")?

because Var%ﬁl = 2a? + a. Therefore by the Assertion 11.1 with § = 5,3, - Eg,%l,
we have

cx (82)] = |ox (82— E82)| = k)2 (2"20) P vardl ke = 3.

Hence - k—2 pt
Cr (XF)| = ®kD? (2"%)" " NVar&} .k = 3,
N 1

because as X2N is a sum of i.i.d. r.v.’s. Apply this and Lemma 11.1 to get: for any
fixed k = 3,4,... and enough large n

)k_z Vargl2

2 k—2
mZNO{(Q‘*‘l) < (k') (2120z) VCH’XIZ,N,

Ck (XF)] = (k)? (2"
since Varg®/2a(a 4+ 1) < 1. Thus r.v. X7 v satisfy the Statulevicius condition

(Sy) withv = 1and A = 213¢, see Appendix. Theorem 11.1 follows from the

Assertion 11.3 with § = X2, 0 = /2Na(a + 1) and A = 213,
Leta < 1.Then

<(2k)'2kZoz < (k) ka < (k)? 21262y g2

p(-em) 28

Hence in this case |Ck ()212\,)| < (kN?*212k-2) Var§12 and |Ck (X%,)| < (k!)?
212Dy rx 12 N-Sorv. X 12  satisfy the Statulevicius condition (S,) with v =
land A = 2!3. Theorem 11.2 follows from the Assertion 11.3 with § =

= /2Na(e + 1) and A = 213,



11 On Tests of Fit Based on Grouped Data 169

Appendix

Let & and &1, &5, ..., beiid. r.v’s with E§ = 0, Varé = 02 > 0, Ci(£) and oy (£)
be respectively, the cumulant and moment of kth order of the r.v. £. The following
two conditions play an essential role in the theory of large deviations (see [35]).
Bernstein’s condition (B,): there exist constants v > 0 and B > 0 such that
lak (6)] < (k)Y FT1B*¥252 forall k =34,....
Statulevicius condition(S,): there exist constants v > 0 and A > 0 such that
ICk(8)] < (k)" A% 52 forall k =3,4,....
Assertion 11.1 ([35]). If £ satisfies condition (B,) then it also satisfies the condi-
tion (Sy) with A = 2B.

Assertion 11.2. Suppose that r.v. £ depending on a parameter A satisfies condition
(S1)G.e., v = 1). Then P {£ > xo} = @(—x) (1 +o(1)) forall x > 0 and x =
0 ((A/o)_l/3) .

Assertion 11.2 is Lemma 2.3 with v = 1 by [35].

Remark. The condition (S,) presented by [35] has the form |Cx(§/0)| < (k!)V*!
A~%=2) then in the Assertions 11.1 and 11.2 one should replace A = ¢/2B by
A=2Bandx = o(AY3) byx = o ((A/U)_l/3), respectively. The slightly
different formulation presented here, of the condition (S,,) is more convenient. For

example, Assertion 11.1 now is easily understood because the following well-known
relation (see, for instance [35, p. 15]).

Assertion 11.3.

k m
_ ar(§) \™
C Zk' -1 my+mo+...+mi—1 Mi4+ms 4+ ... +m 1 1
€)=k (1) (m-+m> - ),Hlml I
where ) _ is summation over all non-negative integer my, ms, . .. , my such that m; +

2my + ...+ kmyp =k
Assertion 11.4 ([31]). For all t: |¢| < 03/n/6B3, with B3 = E |¢]?

] -enl |8l )

E exp
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Chapter 12
Innovation Processes in Logically Constrained
Time Series

Christoph Moller, Svetlozar T. Rachev, Young S. Kim, and Frank J. Fabozzi

Abstract Capturing the relevant aspects of phenomena in an econometric model is
a fine art. When it comes to the innovation process a trade of between a suitable
process and its mathematical implications has to be found.

In many phenomena the likelihood of extreme events plays a crucial role. At
the same time, classical extreme value theory is based on assumptions that cannot
logically be drawn for the phenomenon in question. In this paper, we exemplify the
fitness of tempered stable laws to capture both the probability of extreme events, and
the relevant boundary conditions in a back-coupled system, the German balancing
energy demand.

12.1 Introduction

Classical time series analysis assumes a normal distribution. This assumption is jus-
tified by the central limit theorem (CLT) which states that the infinite sum of random
numbers drawn from any distribution with finite first and second moments con-
verges to the normal distribution. The global financial crisis that began in late 2007,
however, has demonstrated how problematic the assumption of normal innovations
or a Gaussian copular is when it comes to evaluating risk. There is a rich litera-
ture proposing alternative modeling approaches (see, e.g., [21] and the references
within).

In this paper we will analyze a non-financial seasonal autoregressive integrated
moving average (SARIMA) time series whose innovation process is clearly in the
domain of the CLT due to physical boundary conditions. Nonetheless, we will
demonstrate normal innovations to be inadequate in this setting due to the presence
of heavy-tailed distributions in the data. Furthermore, we will address the issue of
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robustness of the Kolmogorow—Smirnow test and the Anderson—Darling test when
testing heavy-tailed distributions.

The remainder of this paper is organized as follows. Section 12.2 reviews clas-
sical linear time series analysis: the non-seasonal autoregressive integrated moving
average (ARIMA) and SARIMA models. Different distribution classes for the corre-
sponding innovation process are discussed in Sect. 12.3 along with distribution tests.
Section 12.4 introduces the balancing energy demand time series and the relevant
boundary conditions. In Sect. 12.5 we describe the SARIMA model used to filter
the innovations that are subsequently analyzed for their distribution in Sect. 12.6.
Section 12.7 concludes and summarizes our results.

12.2 Time Series Models

In econometrics, a linear time series regression can be regarded as a fundamental
building block. We restrict our overview here to the case of discrete time series
models. One widely known example is the autoregressive (AR) model capturing
serial dependence in an analyzed time series. In this model, the realized time series
x; of arandom variable X; is generated from a noise process ¢;, in its standard form
the Gaussian white noise process, and a regression on the past realizations of the
process. One example would be Brownian motion w; in discrete time; however, this
process is not stationary and may grow without limits. Nevertheless, the process is
transformed into a stationary process (w; — w;—_1) by differencing, which in turn
is expanded into the original Brownian motion by summation or integration. So
Brownian motion is a simple example of an integrated stationary AR-process.

If one replaces each past realization in the AR-process by the lagged process, it
is transformed into a possibly infinite regression on past realizations of the noise
process, the so-called moving average (MA) process. Obviously this transformation
is reversible and so any such MA-process can be transformed into a possibly infinite
AR-process. Additionally, Wold’s decomposition theorem states that any stationary
time series may be represented by a possibly infinite MA-process, or equivalently
AR-process. This explains why the combination of the autoregressive moving aver-
age (ARMA)-process has become the standard approach in time series modeling.
The practical advantage over the strict AR- and MA-processes is a parsimonious
usage of parameters. This approach is expanded to an even wider range of non-
stationary time series the ARIMA model, when including the appropriate degree of
differencing to obtain a stationary time series.

Again the consideration of parameter parsimony leads to a further specification,
the SARIMA model. This seasonal ARIMA model is a serial connection of ARIMA
models. Here, one ARIMA model captures a serial dependence structure over cycli-
cal periods (e.g., 1 year), and a further ARIMA model captures the dependence on a
sub-cycle horizon [1]. Using the lag-operator L and the difference-operator V, the
model can be expressed as in (12.1):

dp(L)Pp(L5)VIVPx, = 0,(L)Og(L%)e (12.1)
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Here s is the seasonality, p and P are the orders of the AR-polynomials ¢, @ and ¢
and Q are the orders of MA-polynomials 6 and ®, and d and D are the appropriate
numbers of differencing to obtain stationarity.

Therefore the model is also abbreviated as SARIMA(p,d,q) x (P, D, Q)s.
The crucial difference to the regular ARIMA(p, d, q) model can be best seen by
expanding the multiplicative model into its additive form as in (12.2):

(=1L —poL® — ... = (§s + DL’ — (ps41 — 5P L — . )VIVPy,
=0 4+6,L+6,L%+...4 by + O)LS + (Bs11 + 0,0 LT + . e,
(12.2)

So as an example, a SARIMA(1,0,0) x (1,0,0);, is equivalent to an ARIMA
(13,0, 0) with the constraint ¢13 = —¢d1¢12.

We conclude this section with a brief overview of model identification techniques
that were established by [1]. The first condition a time series needs to fulfill in order
to adapt a linear model is stationarity. This condition is fulfilled if all roots of the
polynomial ¢ (L) lie outside the unit circle. Consequently, the hypothesis of a root
of value one is tested (i.e., the unit root test). Should this hypothesis be rejected, the
time series may be regarded as stationary, otherwise a higher order of differencing
has to be considered. The work of [6] lead to the augmented Dickey—Fuller test
(ADF-test) as an unbiased unit root test.

Once the appropriate degree d of differencing is selected, inspection of the sam-
ple autocorrelation function (SACF) and partial autocorrelation function (SPACF)
will help to select the order of the AR and MA polynomials. The SACF will cut
off at the lag g of a M A(q)-process, while the SPACF will cut off at the lag p of
an AR(p)-process. Unfortunately, the SACF will only gradually tail off for an AR-
process as will the SPACF for a MA-process. Consequently, an A R M A-process will
lead to a tailing off of both SACF and SPACF. Additionally, because it is calculated
on a limited sample, this method is also prone to statistical fluctuations. Nonethe-
less, sudden drops in SACF and SPACF give an indication of appropriate choices
for the parameters p and g of an ARM A-process.

Finally model selection criteria such as the Akaike information criterion (AIC) or
Bayesian information criterion (BIC) should be used as a guide to avoid the pitfalls
of overparameterized modeling.

12.3 Innovations

The main purpose of modeling time series is to identify and understand under-
lying characteristics and then be able to apply this understanding to predict the
future. While a specification of the appropriate parameters of a SARIMA(p, d, q) x
(P, D, Q)s) captures information on the location parameter of a future realiza-
tion, the identification of the distribution of the innovation process ¢; is crucial in
understanding the risk that goes along with anticipating this realization.
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Among all distributions, the class of «-stable distributions stands out as the
distributional class any appropriately scaled sum of independent and identically
distributed (i.i.d.) random variables will converge to. In the context of time series
modeling, this property is of particular interest as the innovations in each time step
are considered to be the sum of many independent events from the same set of
plausible events (i.e., the sum of i.i.d. random variables).

The normal distribution is the most well-known member of the class of a-stable
distributions, and for this distribution the convergence property is known as the CLT.
Relaxing the assumption of existing first and second moments, the CLT is gener-
alized. The limiting distribution is now not generally the normal distribution, but,
instead, a member of the class of «-stable distributions. This is known as the general
central limit theorem (GCLT). In the following we will refer to -stable distributions
as non-Gaussian to distinguish the class from its most well-known member. In gen-
eral, for «-stable distributions there exists no probability density function in closed
form and therefore it is expressed by its characteristic function as given by (12.3):

¢stable(u;a’ O’ /3’ M) = E[eiux] (12.3)
T o
exp (z;m |ou| (1 zﬂ;mgn u) tan > )) a#1
exp (iuu—0|u| (1 + if—(sign u) ln|u|)) ,a =1,
7T

where
1, t>0
signt=4 0, t=0
-1, <0.

In this parameterization, the four parameters (&, 8, o, i) have the following domain
and interpretation:

a: the index of stability or the shape parameter, o € (0, 2)
B: the skewness parameter, f € [—1, +1]

o' the scale parameter, o € (0, +00)

W the location parameter, i € (—o0, +00)

The «-stable distribution is a very rich class of distributions that can capture even
extreme asymmetric and heavy-tailed structural conditions. However, in some prob-
lems the implication of infinite variance, or in the case of « < 1 infinite mean and
variance, is too strong an assumption. At the same time the empirical distribution
might clearly show excess kurtosis or skewness. For these problems the class of tem-
pered stable distributions has been proposed. (See [12,16], and [15]). In an adaption
of the «-stable distribution, the tempered stable distribution has the characteristic
function given by (12.4) with one extra parameter.

pcrs(;a, C Ay, Ao,m) = exp(ium — iuCT (1 —a)(A%1 — 2271 (12.4)
+ CT(—a)(Ay —iw)®* — A% + (A= +iu)® — A%)).
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Here m and C determine the location and scale as do u and ¢ in the a-stable
distribution. However, the skewness is parameterized by A and A_, at the same
time allowing for a faster than «-stable decay in the tails. So a tempered stable
distribution allows for the same flexibility at the center of the distribution as the
«-stable distributions, combined with finite first and second moments. Therefore,
the tempered stable distribution is in the domain of the CLT, though convergence to
the Gaussian limit may be very slow due to the almost «-stable distribution in the
center. So tempered stable distributions are a natural choice whenever the problem
suggests a distribution that is prone to extreme events, but at the same time other
considerations forbid infinite moments; that is, whenever the rate of convergence to
the limiting Gaussian distribution at the infinite sum is too slow to justify a Gaussian
model at the finite time horizon of interest.

The classical tempered stable (CTS) distribution has been introduced under dif-
ferent names in the literature, including the truncated Levy flight by [14], the KoBoL
distribution by [2], and the CGMY distribution by [4].

Having introduced the tempered stable distribution, we conclude this section
with a review of methods to justify the introduction of additional parameters into
a model by specifying a non-Gaussian innovation process. One widely used graph-
ical method is the inspection of the quantile-quantile (QQ)-plot. Figure 12.1 shows
an example of the quantiles of the fitted Gaussian-distribution versus the empiri-
cal quantiles of an observed innovations time series. Ideally, the QQ-plot would be
a straight line of unit slope, and any deviation from this line is an indication of the
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Fig. 12.1 QQ-plot of heavy-tailed data under the Gaussian hypothesis
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Table 12.1 Goodness-off-fit test and their statistic

Test Statistic
KS N1 SUP | Fopp (X) — Fip (x)]
Femp. (X)— Fun.(x)
AD /1 sup ’ o YT Fr 00
o0
CvM n /_OO(FEWIp.(x) - Fth.(x))zdFlhA(x)
00 ( Foup. )= Fu(v) \?
AD? o (m.(’ﬂ(l—m.(n)) dFy.(x)

chosen distribution not adequately describing the innovation time series. One should
keep in mind that because it is based on a finite sample, the empirical quantiles will
never stretch out to arbitrarily small or large quantiles.

Another less subjective method is testing the proposed distribution with a
goodness-of-fit test. All the tests calculate a test statistic (see Table 12.1) to test
the hypothesis of the innovations time series to be a realization of the fitted distri-
bution. These statistics can then be compared to critical values and p-values can be
calculated.

The Kolmogorov—Smirnov (KS) test is known for its critical values to be inde-
pendent of the tested distribution. Like the KS-test, the Anderson—Darling (AD)
test is based on the supremum of the difference of the theoretical and empirical
CDF, however, a weight assigned to each point assigns more weight to the tails of
the distribution. In the case of the «-stable distribution, this feature is obviously of
particular importance.

Both the KS-test and AD-test have to be used with caution when testing a-stable
or tempered stable distributions. We illustrate this drawback in the following exam-
ple. Suppose the underlying distribution is the CTS-(0.3,1,0.5,0.7,0) shown in
Fig. 12.2. There is an almost vertical step in the CDF at the location parameter m.
Because of the dominance of extreme events, the mean of a sample drawn from a
heavy-tailed distribution will only converge slowly to the true mean of zero. Con-
sequently, the steep increase is very likely to be dislocated in the empirical CDF.
In combination with the almost vertical increase at the location parameter, this may
result in large deviances between the empirical and theoretical CDF and a rejection
of the distribution in the KS-test. This argument might even hold true for the AD-test
where the weights are minimal at the location parameter. This behavior is particu-
larly undesirable when analyzing innovation distributions where the true mean of
zero is known and the location parameter is of minor importance. We therefore
suggest a flexibility in adjusting the sample mean before a KS-test or an AD-test.

In contrast to the KS-test and AD-test, the Cramer—von Mises (CvM) test and the
AD?-test are based on the area between the theoretical and empirical CDF (see Table
12.1). The main advantage of these statistics is that they incorporate information
about the total sample, and, in particular, are insensitive to a slight dislocation of the
empirical CDF. The AD?-test introduces a weighting scheme that focuses the test on
the tails of the distribution. One major disadvantage of the CvM-test, AD?-test and
AD-test is that critical values are dependent on the analyzed distribution. One fea-
sible method of obtaining critical values in these cases is to use Monte Carlo-based
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Fig. 12.2 CDF of CTS-(0.3, 1, 0.5, 0.7, 0)

simulation based on the estimated parameter set (see [5]). Therefore, the tests are
not available in common software, and in turn results are less transparent.

12.4 Market Setting and Data

Electricity is a special commodity as it is practically not storable. Consequently, in
electricity grids supply and demand sides have to be in exact equilibrium at all times,
otherwise a blackout will result. This task of active balancing is performed by the
transmission system operator (TSO) who activates bids of up- or down-regulating
energy to maintain equilibrium. In this context, it is important to distinguish between
regulatory energy and balancing energy. Regulatory energy is contracted prior to the
actual balancing action by the TSO to allocate the resources to be able to balance
the grid. Balancing energy is the energy the TSO will settle with balancing respon-
sible entities (BR) who caused a disturbance after the balancing action. The TSO
performs this correction by calling up appropriate regulatory energy bids contracted
prior to the actual balancing.

It is technically not feasible to balance disturbances of arbitrary sizes as the
blackouts in northeast North America in 2003 or the blackout in central Europe
in 2006 demonstrate. So balancing energy demand is constrained around zero;
however, no exact boundary can be specified. In general, balancing energy has to
compensate all unpredictable supply and demand shocks such as a power plant
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outage or a football match going into overtime. Such shocks are unknown in advance
and not predictable over a time horizon of a day or even more. Within the German
electricity market design, balancing energy may however also be used as a substitute
for other energy contracts as long as this position is neither obvious nor exces-
sive (see [3]). This incentive leads to pronounced patterns and serial dependence
(see [19] and [18]).

In this paper we concentrate on the governing innovation distribution of balanc-
ing energy demand in Germany. To uncover this distribution, we therefore have
to filter from the data predictable components. The analytical model used here is
described in [18]. For the German market two factors are identified. These factors
are the gradient of grid load and an arbitrage incentive. Next we restate some of the
results.

The gradient effect is modeled by the out of sample average, which in turn can
be modeled by (12.5) using the load (L(¢)) as a factor.

(VL) = q- (Lq(t) — Lp(t)), ¢ =0.42 (12.5)

This model describes the quarter hourly pattern with a R? of 0.87. The effect is fully
attributable to the discrepancy between the step function of load changes imposed by
the traded contracts on the day-ahead market, and a rather smoothly changing load
in reality. Such effects cannot be observed in most of the other European markets,
because the settlement periods of the day-ahead market and the balancing energy
market are one and the same in these markets (see [9]).

As for the second factor, there exist arbitrage incentives (I(¢), Izec(t)) to sub-
stitute electricity trades on the day-ahead market with strategic positions in the
balancing energy market or avoid technical difficulties in plant operation. This
incentive is driven by the day-ahead prices exceedence of a common price level
and can be modeled as in (12.6). The model captures dependence and saturation
effects, and is compatible with a functional relation that is constant over time as the
parameter estimates and R2-values in Table 12.2 demonstrate.

h(I(t), Lec(t)) = a - ( 2 e~ 1) 4 Liec(t) a € R b,c € Ry (12.6)

1+b-e

Table 12.2 Parameters and R? fitting to out of sample data. Source [18]

Parameters R? factor model
Year a[MWh] b[E] c[€7'] I(t)only I(t)and I,..(t)
2003  940.045 1.053  0.035 0.6948 0.7252
2004 901.082 1.113  0.039 0.4448 0.6170
2005 918.633  1.089  0.039 0.6069 0.7518
2006 902.485 1.072  0.038 0.8424 0.8499
2007  928.798 1.081  0.037 0.7571 0.7998

2008  884.110 1.068  0.043 0.6530 0.7025
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The sample variance is reduced by 12 and 19% in the case of the gradient effect
and the arbitrage incentive, respectively. This clearly demonstrates the existence of
non-random predictable components in the balancing energy demand data.

The raw data are obtained from the publications of balancing energy demand
the four German TSOs from January 2003 to December 2008. Namely the four
sources are RWE [22], E.on [8], EnBW [7], and Vattenfall [24]. These data from
four sub-zones are combined to obtain one hypothetical German zone to account
for interzonal balancing effects and provide a better correspondence to the single
German day-ahead market EEX.

12.5 SARIMA Model of Balancing Energy Demand

Here we focus on the analysis of the residual time series after subtracting the effects
described by the analytical model given by (12.5) and (12.6). Before trying to adapt
a linear time series model to the residuals, the data need to be checked for station-
arity. We apply an augmented Dickey—Fuller unit root test. The null hypothesis of
a unit root is rejected at a significance level below o = 0.001 even when including
the first 168 lags for the regression.

This finding is supported also by a consideration of the physical boundary con-
ditions of the underlying data. Balancing energy demand is fulfilled by the grid
operator to ensure grid balance. This energy has to be delivered physically by power
stations, and so the installed capacity imposes a hard boundary. This boundary can,
however, never be reached, as the response time and response capacity of power sta-
tions impose an even tighter boundary. Due to their design, powers stations cannot
run on an arbitrary fraction of their designed capacity, but instead have to be oper-
ated within a certain bandwidth. Additionally, a complex system such as a power
station has a considerable amount of inertia, and cannot instantaneously adapt to
changes in operation. When looking at the total generation stock, although these
facts do not translate into a hard boundary, the true limits will depend on the exact
condition and history of all individual facilities connected to the grid. Nonetheless a
limit to fluctuations the grid operator can manage always exists. So it is physically
impossible for the balancing energy demand to grow to very large positive values
or fall to very small negative values, but balancing energy will always be within a
bandwidth around zero. Mathematically, this argument relates to a stationary time
series, and the absence of unit roots. We can therefore model the data without the
need of further differencing.

As a first step, an inspection of the SACF and SPACF of the residuals in Fig. 12.3
shows the presence of SARIMA effects in the data. The autocorrelation decays off
with increasing lag. Additionally, this decay is disturbed at multiples of 24, indi-
cating a seasonality of 24. This picture is supported by the partial autocorrelation
function displaying a drop at lag one and 24, together with a decaying negative
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Fig. 12.3 Sample autocorrelation and partial autocorrelation

Table 12.3 Parameter estimates of SARIMA model

SARIMA
(1,0,0) X (1,0, 1)24 (3,0,0) X (1,0, 1)24
Gaussian t(v) Gaussian t(v)
ary 0.8185 0.8238 0.7974 0.8036
ar; - - 0.0084 0.0079
ary 0.9572 0.9571 0.9419 0.9427
mayy —0.8502 —0.8529 —0.8291 —0.8328
o 341.9658 341.9410 341.7744 342.0834
v - 9.1080 - 9.1396
AIC 763,170 762,080 763,060 761,970
BIC 763,200 762,110 763,090 762,000

partial autocorrelation at lags following multiples of 24. Moreover, the SPACF indi-
cates another step at lag three. We therefore choose SARIMA(1,0,0) x (1,0, 1)24
and SARIMA(3,0,0) x (1,0, 1)24 as candidates for the model. Additional to the
classical model with Gaussian innovations we include models with ¢-distributed
innovations as representatives of heavy-tailed innovations in the analysis. This
approach is suggested by [25] as a compromise between a heavy-tailed innova-
tion distribution and robust parameter estimates for the SARIMA-model. Table 12.3
holds the AIC and BIC values of different specifications, including both Gaussian
and 7-distributed innovations. We choose the SARIMA(1, 0, 0) x (1,0, 1)24 model
with ¢-distributed innovations for two reasons. First, the AIC and BIC values indi-
cate a preference of ¢-distributed innovations over the Gaussian case. Second, the
ars coefficient is small and, as we will discuss below, coefficients at low lags are
of minor practical relevance. Note that the 7-distributed innovations demonstrate the
necessity of a heavy-tailed noise term in the model. This will be further investigated
in Sect. 12.6.

We conclude this section with an analysis of the consistency of the model over
time and a test of its forecasts. Table 12.4 reports the parameter estimates of the
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Table 12.4 Parameter estimates of SARIMA model

Parameter a ayy by o v
Total 0.8238 09571 —0.8529 341.9410 9.1080
2003 0.8522 0.9307 —0.7973 341.9410 9.4983
2004 0.7860 0.9421 —0.8184 328.8776 12.3724
2005 0.7810 09359 —0.8248 325.6431 10.7623
2006 0.8092 0.9455 —0.8397 336.8019 9.0538
2007 0.8067 09586 —0.8620 329.9785 10.1529
2008 0.7861 09515 —0.8541 341.9410 9.2676

model based on yearly sub-samples. The parameter estimates are very consistent
with the overall model. We therefore decided to test the forecasts of the overall
model rather than the individual yearly models.

The information on balancing energy demand is not continuously revealed to the
market, but rather published only once a month, including the data for the preceding
month. So the data for May will be available by July. We therefore do not test the
one-time step forecast, as this has no practical implication in this market. Instead,
we test a forecast adapted to the information revealed to the market. As aresult, fore-
casting is performed once a month based on the information lagged 1 month (i.e.,
the forecast horizon is 720-1,440 lags). Because one of the TSOs also publishes the
balancing demand data in its zone with a time delay of only 3 days, we also test
the implications of the model on this time horizon (i.e., a forecast horizon of 72-96
lags). In both cases that we test, the sample variance is reduced by subtracting the
conditional expectation. When using the monthly forecast, the variance is reduced
by 3.69%; applying a 3-day forecast horizon results in a 11.22% reduction.

This additional variance reduction as compared to the analytical model in
Sect. 12.4 can be decomposed into two components. For the first component there
exist comparatively short-lived patterns in the data. These patterns can be under-
stood as a linear correction term for the analytical model. The second component
captures a non-zero conditional mean of the time series. Neither the gradient effect
nor the arbitrage incentive described in Sect. 12.4 can explain a non-zero expectation
value of the balancing energy demand over extended periods of time. Both effects
will average to zero over a few cycles of their respective seasonality. However when
looking at the average forecast of the SARIMA-model in Table 12.5, it is evident
the SARIMA forecast does not average to zero over a few cycles. Furthermore, it
is argued in [17] that this non-zero mean over extended periods of time is sufficient
to influence the electricity price on the day-ahead market. Other studies of market
power abuse in Germany identify the same periods as periods of abuse, which show
highly negative mean SARIMA forecasts (i.e., artificial demand) in their analysis
(see [10,11,23]).
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Table 12.5 Average prediction of SARIMA model
Average prediction

Horizon 2003 2004 2005 2006 2007 2008

One month  —198.9537  —179.9403 —110.6992  21.8594 —118.4479  —52.1579
Three days  —372.3728  —307.2270 —214.6126  18.6222 —217.6485 —190.4806

12.6 Innovation Distribution

The parameter estimation of the SARIMA-model used in Sect. 12.5 is based on a ¢-
distributed innovation process. Models with Gaussian innovations were disregarded
based on AIC and BIC values reported in Sect. 12.5. In this section, we will further
investigate the heavy-tailed innovation process.

The innovations process is of particular importance in the balancing energy mar-
ket as it governs the risk involved with balancing the grid. In general, TSOs have to
allocate sufficient regulatory power to be able to maintain grid operation and avoid a
blackout. The capacity that is considered sufficient is usually defined by a threshold
probability for a blackout (i.e., the probability of fluctuations exceeding the allo-
cated capacity). So the more precise the quantiles of the innovations distributions
are known, the more efficiently resources may be allocated.

The first step of the investigation is the QQ-plot of the innovations time series
and the fitted -distribution in Fig. 12.4. From this figure it can be seen that the
t-distribution does not adequately capture the risk in the tails of the empirical
distribution because the QQ-plot deviates from the diagonal.

Due to the conceptual advantage of modeling the data with a distribution in the
proximity of the GCLT as discussed in Sect. 12.3, we test both the «-stable and the
CTS-distribution as a more adequate model for the innovations time series. Both
distributions are estimated by the Fourier inversion formula and their characteristic
functions given by (12.3) and (12.4). This inversion is, in turn, numerically esti-
mated by the fast Fourier transform (FFT) method. A more detailed description of
the method is given in [20] and [13]. Table 12.6 shows the estimated parameter sets.
As can be seen in Fig. 12.5, the heavy-tailed distribution captures the likelihood of
extreme events more accurately than the ¢-distribution.

In the next step, all three distributions are compared using the goodness-of-fit
tests mentioned in Sect. 12.3. The results are summarized in Table 12.7.

The p-values of the KS-test clearly indicate that the CTS-distribution describes
the innovations best, as its p-value is 49 and 9 orders of magnitude greater than
the p-value of f-distribution and «-stable distribution, respectively. However the
p-value of the CTS-distribution is still low. As discussed in Sect. 12.3, the KS-test
is responsive to small fluctuations in the location parameter, while such fluctuations
are to be expected with heavy-tailed distributions. Also, the SARIMA model implies
a location parameter of zero for the innovation process, so we do not need to focus
on the location parameter. We therefore correct the mean of the innovations time
series for such fluctuations within the 95% confidence bounds. The corresponding
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Fig. 12.4 QQ-plot ¢-distribution

Table 12.6 Estimated parameters of heavy-tailed distributions

Distribution Parameters

«-stable « o p ®
1.9107 0.0048 0.6711 0-fixed

CTS o C Ag A m
0.9122 1.4856 1.5168  O-fixed

[ —
Ie—a)(A242072)

statistics are identified by an asterisk (*). Again the CTS-distribution provides the
best description of the data. Furthermore, the CTS-distribution is acceptable at a 5%
significance level. The other statistics reported provide further support for selecting
the CTS-distribution over both the ¢-distribution and the «-stable distribution.

12.7 Conclusion

The wide application of linear time series models in finance has made the short-
comings of inadequate innovation processes and correlation structures evident.
Tempered stable distributions have been proposed and tested on financial time series
data to overcome these problems. The tempered stable distribution is an expan-
sion of the a-stable distribution. It combines heavy-tailed innovations over multiple
timescales with finite higher moments. These properties make tempered stable
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Table 12.7 Goodness-of-fit statistics and p-values
Distribution
t a-stable CTS
Test Statistic p-value Statistic p-value Statistic p-value
KS 0.0331 1.37-10—% 0.0156 1.75-10~1 0.0082 0.0016
AD 0.0883 - 0.0333 - 0.0170 -
KS* 0.0316 5731074 0.0134 1.40-107% 0.0059 0.0538
AD* 0.0854 - 0.0316 - 0.0126 -
CvM* 23.4809 - 2.2939 - 0.5144 -
AD?x 625.3888 - 685.0225 - 611.5575 -

distributions an excellent choice for modeling phenomena dependant on extreme
events, which are at the same time bounded by other considerations.

In this paper, we apply the classical tempered stable model to German balanc-
ing energy demand data and demonstrate its fitness. The CTS-distribution describes
the risk of unpredictable events in the electricity grid, while at the same time cap-
turing physical boundary conditions in the model. In the balancing energy market,
these advantages can be used to allocate the resource capacity more efficiently, and
thereby reduce grid tariffs. Additionally, the SARMIA model is able to separate
predictable balancing energy demand from unpredictable shocks. In general, the
predictable fraction of balancing energy demand can be satisfied by a wider and
technically less demanding range of providers. The German market design includes



12 Innovation Processes in Logically Constrained Time Series 187

the option to trade this predictable fraction in strategic positions on the balancing
market, while the reserve capacity for the unpredictable shocks is traded on the
market for regulatory energy. A further development of strategic positions in the bal-
ancing energy market could therefore increase the competitiveness of the capacity
reserve market. In view of the planned increase of highly fluctuating regenerative
power in the German system, this advantage will become even more pronounced in
the future.
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Chapter 13
Laws of Large Numbers and Nearest Neighbor
Distances

Mathew D. Penrose and J.E. Yukich

Abstract We consider the sum of power weighted nearest neighbor distances in a
sample of size n from a multivariate density f of possibly unbounded support. We
give various criteria guaranteeing that this sum satisfies a law of large numbers for
large n, correcting some inaccuracies in the literature on the way. Motivation comes
partly from the problem of consistent estimation of certain entropies of f.

13.1 Introduction

Nearest-neighbor statistics on multidimensional data are of long-standing and con-
tinuing interest, because of their uses, e.g., in density estimation and goodness-of
fit testing [3, 11,21], and entropy estimation [2, 4,9, 10]. They form a multivariate
analog to the one-dimensional spacings statistics in which the work of S.R. Jam-
malamadaka, the dedicatee of this paper, has featured prominently. For example,
[16] uses nearest neighbor balls to generalize the maximum spacings method to
high dimensions and to establish consistency in estimation questions.

In the present note we revisit, extend and correct some of the laws of large num-
bers concerned with sums of power-weighted nearest-neighbor distances that have
appeared in recent papers, notably [10, 15, 19].

Fix d € Nand j € N. Given a finite X C R4, and given a point x € X, let
card(X) denote the number of elements of X', and let D(x, X) := D;(x, X) denote
the Euclidean distance from x to its jth nearest neighbor in the point set X' \ {x},
if card(X) > j; set D(x, X) := 0if card(X) < j. Let f be a probability density
function on R¥, and let (Xi)ien be a sequence of independent random d -vectors
with common density f. Forn € N, let X, := {X1,..., X,}. Let € R and set
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Snai= Y D0, %)% =) (DX, X))

XEXy i=1

Certain transformations of the S, o have been proposed [9, 10] as estimators for
certain entropies of the density f which are defined in terms of the integrals

= [ rordy (>0,

For p > 0 with p # 1, the Tsallis p-entropy (or Havrda and Charvit p-entropy
[7]) of the density f is defined by H,(f) := (1 —1,(f))/(1 — p), while the Rényi
entropy [17] of f is defined by H(f) :=log I,(f)/(1 — p).

Rényi and Tsallis entropies figure in various scientific disciplines, being used in
dimension estimation and the study of nonlinear Fokker—Planck equations, frac-
tal random walks, parameter estimation in semi-parametric modeling, and data
compression (see [4] and [10] for further details and references).

A problem of interest is to estimate the Rényi and Tsallis entropies, or equiv-
alently, the integrals I,(f), given only the sample {X;}7_, and their pairwise

distances. Let wg = 7%/2/I'(1 + d/2) denote the volume of the unit radius
Euclidean ball in d dimensions, and set y(d, j) := a);“/ d (W) This note

provides sufficient conditions on the density f establishing that y(d, j) ™ 'n"1S, o
converges t0 I1_q/4(f) in L, or in L2, In other words, since L' convergence
implies convergence of means, we provide sufficient conditions on f guaranteeing
that y(d, j)"'n"18, 4 is an asymptotically unbiased and consistent estimator of

Ii—gra(f).

13.2 Results

Two of our results can be stated without further ado.

Theorem 13.1. Let o > 0. Suppose the support of f is a finite union of convex
bounded sets with nonempty interior, and f is bounded away from zero and infinity
on its support. Then as n — oo we have L? and almost sure convergence

als mald (F(j +a/d)
“ Q)

Theorem 13.2. Letq = 1 orq = 2. Let o € (—d /g, 0) and suppose f is bounded.
Then (13.1) holds with L4 convergence.

)Il—a/d(f)- (13.1)

For the interesting case when o > 0 and f has unbounded support, our results
require further notation. Let | - | denote the Euclidean norm on R<. For r > 0, define
the integral
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M) =EX] = [l o,

and define the critical moment r.( f) € [0, co], by

re(f) :=sup{r >0: M,(f) < oo}.

If r < s and Ms(f) < oo, then M,(f) < oo. Hence M, (f) < oo forr < r.(f)
and M, (f) = oo forr > r.(f).

For k € N, let A; denote the annular shell centered around the origin of R4
with inner radius 2% and outer radius 2"“, and let Ay be the ball centered at the
origin with radius 2. For Borel measurable A C R¥, set F(A) := P[X, € A] =
J4 f(x)dx.

We can now state the rest of our results.

Theorem 13.3. Letq = 1 0rq = 2. Leta € (0,d/q). Suppose I1_yq(f) < 0o,
andre(f) > qad/(d — qa). Then (13.1) holds with L4 convergence.

We shall deduce from Theorem 13.3, that when f(x) decays as a power of |x|,
the condition /;_q/4(f) < oo is sufficient for L' convergence:

Corollary 13.1. Suppose there exists B > d such that f(x) = O(|x|™#) as |x| —
00, i.e., such that for some finite positive C we have

CUxI™ < f(x) <C|x|™, VxeR?, |x|=C. (13.2)

Suppose also that I1_g/q(f) < oo for some a € (0,d). Then (13.1) holds with L'
convergence.

Our final result shows that in general, the condition /1_4/4(f) < oo is not
sufficient alone for L! convergence, or even for convergence of expectations. It can
also be viewed as a partial converse to Theorem 13.3 showing, under the additional
regularity condition (13.3), that when ¢ = 1 the condition r.( ) > qad /(d — qa)
is close to being sharp.

Theorem 13.4. Let0 < o < d. Then (i) If rc(f) < ad/(d — ), and also for some
ko € N we have

. F(Ax) F(Ax)
0< inf ——— < sup ——— < o0, (13.3)
k>ko F(Ag-1) ~ k>ko F(Ak-1)

then lim sup,,_, oo E[n71S, o] = 00;

(ii) For 0 < r < ad/(d — a) there exists a bounded continuous density function
f on R? satisfying (13.3), such that Ii_g/a(f) < oo, but with re(f) = r so that
limsup,,_, o E [n71 Sy o] = 00 by part (i).

The value of the limit in (13.1) was already known (see Lemma 13.1). The con-
tribution of the present paper is concerned with the conditions under which the
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convergence (13.1) holds; in what follows we compare our conditions with the exist-
ing ones in the literature and also comment on related limit results. For conditions
under which n=1/2 (Sn,a —E Sy o) is asymptotically Gaussian, we refer to [1,12,14].

Remark. (i) Theorem 13.1. The condition in Theorem 13.1 is a slight relaxation of
condition C1 of the L? convergence results in [15] or [19], which assume a poly-
hedral support set. When the support of f is the unit cube, Theorem 2.2 of [8]
gives an alternative proof of almost sure convergence in (13.1) (we remark that
Theorem 2.2 of [8] contains an extraneous [E in the left-hand side). Theorem 8.3
of [20] uses subadditivity to obtain (13.1) when the support of f is the unit cube
and when o = 1, but it does not identify the limiting constant. The convergence
of means implied by Theorem 13.1 was previously obtained, under some extra
differentiability conditions on f, in [5].

(ii) Theorem 13.2. The L' convergence of Theorem 13.2 improves upon Theorem
3.1 of [10], which establishes mean convergence; the L? convergence of Theorem
13.2 is contained in Theorem 3.2 of [10] and we include this for completeness.

(iii) Theorem 13.3. The condition in Theorem 13.3 corrects the condition of the
corresponding result given [15], where for L' convergence it is stated that we need
re(f) > d/(d — a); in fact we need instead the condition r.(f) > ad/(d — ).
In the proof of Theorem 13.3 below, we shall indicate the errors in the proof in
[15] giving rise to this discrepancy. This correction also applies to condition C2 in
Theorem 2 of [19], the proof of which relies on the result stated in [15].

(iv) Theorem 13.4. The condition (13.3) holds, e.g., if f(x) is a regularly varying
function of |x|. Given (13.3) and given /;_q/q < o0, Theorem 13.4 shows that
the condition r.(f) > ad/(d — &) is necessary for L' convergence of n™1S, 4,
while Theorem 13.3 says that r.(f) > ad/(d — «) is sufficient. It would be of
interest to try to find more refined necessary and sufficient conditions when r.( ) =
ad/(d —a).

(v) General ¢. For ¢ : Rt — R* put S, 4 = ZXEX}'! ¢4 D(x, X,)). If
¢ has polynomial growth of order «, that is if there is a constant & € (0, co) such
that ¢(x) < C(1 4+ x%) for all x € RT, then straightforward modifications of the
proofs show that under the conditions of Theorem 13.1 or Theorem 13.3 we have
the corresponding L4 convergence

7 Sup > [ EDDO.Psco) (i

where for all z > 0, P, is a homogeneous Poisson point process in R? having
constant intensity t, and D(0, P;) is the distance between the origin of R4 and its
Jj th nearest neighbor in P;.

(vi) Minimal spanning trees. Given a finite ¥ C R? and ¢ : RT — R, let

Ly(X):= D (e,

eeMST(X)
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where MST(X) denotes the edges in the graph of the minimal spanning tree on X’
Thus Lg(X) is the sum of the ¢-weighted edge lengths in the minimal spanning tree
on X.Letg = 1 or 2. If ¢ has polynomial growth of order «, with @ € (0,d/q), if
Ii—qja(f) <oo,andif re(f) > qad/(d — qo) then, as may be seen by following
the proof of Theorem 13.3, the proof of Theorem 2.3(iii) of [15] in fact shows that
as n — oo we have

1
Loty =5 [ E| X aled | st

R4
e€MST(0,P r(x))

where the convergence is in L4, and where MST (0, P f(x)) denotes the edges in the
minimal spanning tree graph on 0 U P r(y) incident to 0, the origin of R?. When
q = 2, this is new whereas for¢ = 1 and @ € (0, 1), this improves upon Theorem
2.3(iii) of [15], which requires r.( f) > max(ad/(d — @), d/(d — )).

(vii) Non-existence of density. If the {X;}"_, fail to have a density, then nor-
malization of S, , may involve exotic functions of n, including log periodic nor-
malizations, as is the case when the {X;}”_, have a Cantor distribution on [0, 1];
see [18].

(viii) Comparison with [10]. The convergence of expectations corresponding
to (13.1) is given as the main conclusion in Theorem 3.1 of [10]. In the case
1 —a/d < 1 of that result, it is claimed that this convergence of expectations
holds without any extra conditions besides finiteness of I;_q /4. Theorem 13.4 here
disproves this assertion; the argument in [10] requires that convergence in distribu-
tion implies convergence of rth moments, which is not in general true. On the other
hand, Corollary 13.1 shows that if we assume f(x) decays as some power of |x|
then finiteness of /;_y/4 is indeed a sufficient condition for convergence in L', and
hence also convergence of expectations.

13.3 Proofs

This section provides the proofs of the results stated in the preceding section. We
denote by ¢, C,C’, and C” various strictly positive finite constants whose values
may change from line to line. The proofs of Theorems 13.1, 13.2 and 13.3 use the
following result.

Lemma 13.1. Let g € {1,2} and o € R. Suppose for some p > q that E[(n'/¢
D (X1, X,))*P] is a bounded function of n. Then (13.1) holds with L9 convergence.

Proof. Since D is a stabilizing functional on homogeneous Poisson point processes
[15], we can apply Theorem 2.2 of [15] or Theorem 2.1 of [15] to get LY conver-
gence of n~1S, 4 to a limit which is expressed as an integrated expectation in [15]
(see (2.15) of [15]). It was shown in [19] that this limit is equal to the right hand
side of (13.1) (and this is also consistent with the limiting constant in [5]). O
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Proof of Theorem 13.1. Recall that we assume the support of f, namely supp
(f) == {x € R? : f(x) > 0}, is a finite union of bounded convex sets
with nonempty interior, here denoted Bji,..., By,. Set A := sup{|x — y| : x €
supp(f), y € supp(f)}, the diameter of the support of f. By assumption, A < oo.
Also we assert that there is a constant ¢ > 0 such that for r € (0, 4],

F(Br(x)) > cr?, Vx e supp(f). (13.1)

To see this, take §; > 0 such that for 1 < i < m there is a ball B;” of radius §;
contained in B;. There is a constant 55 > 0 such that for 1 <i < m, if x € B;, and
r < 81, then the intersection of the ball of radius r centered at x with the convex
hull of the union of B;” and x has volume at least 52rd . This region is contained
in B; and (13.1) follows for r € (0, §1]. But then (with a different choice of ¢)
(13.1) follows for r < A. Hence, for 0 < ¢t < An'/4 and with B(x;r) denoting the
Euclidean ball of radius r centered at x,

Pn"iD(X,,X,) >t] < sup Pleard(X,—; N B(x;n" V1)) < j]

xesupp(f)
i n—1
< _ (cn_ltd)i(l _ Cn—ltd)n—l—l'
(%)
j—1
<C Ztid exp(—cn_ltd(n —1-1))
i=0

< C'(1 + 1Y) exp(—ct?) < C" exp(—(c/2)t%).

Moreover this probability is clearly zero for t > An'/?. Hence, for & > 0 and
p>2,

o0
B[ D)) = [P DG ) > Pl
0
o0
<C [ exp(—(c/2)u’@P)yay
0

which is finite and does not depend on n. Therefore we can apply Lemma 13.1 to
get the L2 convergence (13.1).

For almost sure convergence, we apply Theorem 2.2 of [13], where here the test
function considered in that result (and denoted f there, not to be confused with
the notation f as used here) is the identity function. It is well known (see [3], or
Lemma 8.4 of [20]) that there is a constant C := C(d) such that for any finite
X C R4, any point x € X is the jth nearest neighbor of at most C other points
of X. Therefore adding one point to a set X within the bounded region supp( /)
changes the sum of the power-weighted j th nearest neighbor distances by at most a
constant. Therefore (2.9) of [13] holds here (with § = 1 and p’ = 5 say), and the
almost sure convergence follows by Theorem 2.2 of [13]. O
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Proof of Theorem 13.2. The proof depends on the following lemma. Recall that
(X;)i>1 are ii.d. with density f. Given X;, let V, denote the volume of the
d -dimensional ball centered at n'/4 X 1 whose radius equals the distance to the jth
nearest point in n'/¢ (X, \ X;), where for r > 0 and X C R¢ we write r X for
{rx:x € X}.Forall x € R4, foralln =2.3,...and forall v € (0, 00) let

Fux(v) = PV, <v|X1 =x]. (13.2)

Lemma 13.2. If f is bounded and § € (0, 1), then
o0
supE Vn_5 = sup/ [ v_‘ngn,x(v)f(x)dx < 00.
n n JrRA Jo

Proof. Since [;° v PdF(v) = p [ v P~ F(v)dv for any p € (0,1) whenever
both integrals exist (see, e.g., Lemma 1 on p. 150 of [6]), we have for all x € R4

o0 o0
/ v—ﬁdF,,,x(v)zsf VIR, c(dy
0 0
1 oo
f/ v_s_an,x(v)dv—l—S/ v 1y
0 1
1
:/ VIR, c(dy + 1.
0

With B, (x) denoting the ball of volume v around x, for all v € (0, 1) we have

Fax(v) = P[Va vIX1 = x] = 1 = Pleard(n'/* %, 1 B,(n"/x) < j]
1 — Plcard(n'/? X, N B,(n'/9x)) = 0]

n—1
1_(1_ / f(z)dz) . (13.3)
B,/n(x)

Since f is assumed bounded we have

A

Fnx(v) = 1—exp ((n — 1) log(l — || f loov/7)) .

When n is large enough, for all v € (0, 1) we have (n — 1) log(1 — || f|leov/n) >
—2| f looV, and so for all x € R?

Fox(v) = 1T =exp(=2[ flloov) = 2/ f lloov.

Hence for all n large enough and all x we have fol VvIITIE,  Mdv < 2| flleo

fol v8-1yq v, demonstrating Lemma 13.2.
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Now to prove Theorem 13.2, we choose p > ¢ such that —1 < ap/d < 0 and

invoke Lemma 13.2 to conclude sup,, E [V;;"? / d] < 00. We now apply Lemma 13.1
to complete the proof of L? convergence. O

The proof of Theorem 13.3 uses the following lemma. Recall from Sect. 13.2 the
definition of the regions A,k > 0.

Lemmal3.3. Let 0 < s < d. If re(f) > sd/(d — s), then Y 5o, 2k
(F(Ap))@=9/d < .

Proof. We modify some of the arguments on p. 85 of [20]. For all ¢ > 0, by Holder’s
inequality we have

Z 2ks (F[Ak])(d—s)/d — Z 2—£ks (F[Ak])(d—s)/d2(l+8)ks
k k

s/d
< (Z(z—ekS)d/S) (Z F[Ak](z(l-‘re)kS)d/(d—s))
k k

(d—s)/d

(d—s)/d
< C(e.s) (Z fA |x|“+6>~‘d/<d—~‘>f<x>dx)
k k

which, for ¢ small enough, is finite by hypothesis. O

Proof of Theorem 13.3. We follow the proof in [15], but correct it in some places
and give more details in others. We aim to use Lemma 13.1. Since we assume 0 <
a < d/q,we can take p > g with ap < d. Clearly

E[(n"4D(X;, X,))*?] = n®P/471E [Z D(X,-,Xn)"‘p:| = n®P/TIE [LP (X)),
i=1

(13.4)

where for any finite point set X C R?, and any b > 0, we write L?(X)
for D v D(x, X)? (and set L?(¥) := 0). Note that for some finite constant
C = C(d, j) the functional X — L?(X) satisfies the simple subadditivity relation

LP(xuY) < LP(x) + L) + Cct® (13.5)

for all # > 0 and all finite X and ) contained in [0, t]d (cf. (2.2) of [20]).
Asin (7.21) of [20] or (2.21) of [15] we have that

oo

L% (X,) < <§ LP(X N Ak)) +C(p) max X, (136)
<i<n
k=0 -

In the last sentence of the proof of Theorem 2.4 of [15] it is asserted that the last
term in (13.6) is not needed, based on a further assertion that one can take C = 0
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in (13.5) here, but these assertions are incorrect. For example, if card())) < j then
LP(Y) = 0 but L2(X U Y) could be strictly greater than L?(X). Similarly, if
card(X;,, N Ax) < j then the term in (13.6) from that k is zero but the corresponding
contribution to the left side of (13.6) is non-zero.

Combining (13.6) with (13.4) yields

E[(n'/? D(X1, X,)*?] < n@r=DI9E [Z L7 (X, 0 Ak)}
k
+ C(p)E [n©@P=D/4 max | X;|*P]. (13.7)
1

By Jensen’s inequality and the growth bounds L*P(X) < C(diamX)*?
(card(X))@—eP)/d (see Lemma 3.3 of [20]), we can bound the first term in the
right hand side of (13.7) by

szkap(F[Ak])(d—Otp)/d_ (13.8)
k

Recall that we are assuming 0 < & < d/q and also r.(f) > gda/(d —qa) (the last
assumption did not feature in [15], but in fact we do need it). Let p > g be chosen
sothatre(f) > dap/(d —ap) as well as ap < d. Setting s = ap in Lemma 13.3,
we get that the expression (13.8) is finite. Thus the first term in the right hand side
of (13.7) is bounded by a constant independent of n.

The second term in the right hand side of (13.7) is bounded by

1
C(p) ([ g |: max |X;|*? > tn(d_ap)/di| dt
0

1<i<n

o
+/ P |:max | X; %P ztn(d_“p)/di| dt)
1

1<i<n

o0
<C(p) (1 +n/1 P|X, [#Pd/(@=ap) > ,d/(d—ap)n]d,).

By Markov’s inequality together with the assumption r.(f) > dap/(d — ap), this
last integral is bounded by a constant independent of n.

Therefore the expression (13.7) is bounded independently of n, so we can apply
Lemma 13.1 to get the LY convergence in (13.1). O

Proof of Corollary 13.1. Suppose for some 8 > d that f(x) = O(|x|7#) as |x| —
oo. Then it is easily verified that given a € (0, d), the condition /;_q/q(f) < 00
implies that —(1 — «/d) + d < 0 and hence B > d?(d — a)~'. Moreover,
it is also easily checked that r.(f) = B — d so thatif 8 > d?(d — )~ then
re(f) > da/(d —a).

Therefore, if I1_4/4(f) < 0o we can apply the case ¢ = 1 of Theorem 13.3 to
get (13.1) with L! convergence. O
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The proof of Theorem 13.3 shows that

supE [(n"? D(X1, X,))?] < 0. (13.9)

if ¢ > Ois such that ed /(d — &) < rc(f). The proof of Theorem 13.4, given below,
shows that the condition ed /(d — €) < r.(f) cannot be dropped in general.

Proof of Theorem 13.4. Let 0 < @ < d. Suppose that r.(f) < ad/(d — «), and
(13.3) holds for some k¢ € N. Choose r,s such that r.(f) <r < s < ad/(d —
@). Then M, (f) = o0, 50 ) 2"k F(Ay) = oo and therefore there is an infinite
subsequence /C of N such that

2k F(A) =1, kek. (13.10)

Indeed, if no such KC existed, then for all but finitely many k we would have
27"k F(Ag) < 209k which is summable in k.

Given k € N, and set n(k) = [(F(Ax))~'], the smallest integer not less than
(F(Ag))™!. Let E be the event that X; € Ay but X; ¢ Ax_; U Ay U Apy; for
2 <i < n(k). Then by the condition (13.3), there is a strictly positive constant c,
independent of k, such that for k > ko we have

P[Ex] = F(Ax)(1 = F(Ag—1 U A U A 11))"®71 > cF(A4p).

If Ej occurs then D(X1, X)) > 2k=1 5o for n = n(k) we have (for a different
constant ¢) that

E[n~'Spa] = E[(nY9D(X1, X,))%] = n¥9E[D(X1, X,)*1E, ]

> en®4F(Ar)2 = c(F(Ap))' ™92k,
By (13.10), for k € I this lower bound is at least a constant times 2k(a—s(d—a)/d)
and therefore tends to infinity as k — oo through the sequence /C, concluding the
proof of part ().

For part (ii), for each k > 2 choose, in an arbitrary way, a unit radius ball By
that is contained in Ag. Given r € (0, ad /(d —)), consider the density function f
with f(x) = C27"* for x € B,k > 2, and with f(x) = 0 forx € R? \ U By;
here the normalizing constant C is chosen to make f a probability density function.
This gives F(Ax) = Cwgy27 "% for each k > 2; it is easy to see that this f has
r¢(f) = r, and that (13.3) holds with kg = 3. Also, forany p > O we have I,(f) =
®gCP Y 1on 277Pk which is finite, so in particular /;_, /a < oo. This choice of f is
bounded but not continuous, but can easily be modified to a continuous density with
the same properties, e.g., by modifying f in an annulus near the boundary of each
ball By so as to make it continuous, and then adjusting the normalizing constant C
accordingly. O
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Chapter 14

Nonparametric and Probabilistic Classification
Using NN-balls with Environmental

and Remote Sensing Applications

Bo Ranneby and Jun Yu

Abstract National and international policies today require environmental follow-
up systems that detect, in a quality assured way, changes over time in land use and
landscape indicators. Questions related to environmental health and spatial patterns
call for new statistical tools. We present in this paper some new developments on the
classification of land use by using multispectral and multitemporal satellite images,
based on techniques of nearest neighbour balls. The probabilistic classifiers intro-
duced are useful for measuring uncertainty at pixel level and obtaining reliable area
estimates locally. Also some theoretical considerations for the reference sample plot
method (today named k-NN method in natural resource applications) are presented.

14.1 Introduction

Environmental pressures on ecosystems and the continuous loss of suitable habi-
tats for different species jeopardize the ambitions of a sustainable development.
Environmental work needs to be enforced at a global, national, regional and local
level.

Spatial patterns at different scales are important characteristics of environmental
quality. Spatial elements that indicate high environmental quality are usually quite
rare and important changes are sometimes very subtle. Existing monitoring pro-
grams linked to landscape surveillance do not permit evaluation of data at a local
level or of smaller regions. The landscape perspective requires wall-to-wall analysis
that are scale-independent, i.e., the results can be used at different levels and the
accuracy of the measurements can be clearly defined.

Remote sensing of satellite images offers great potential to assess wall-to-wall
changes in the health of ecosystems and identify risks. So far, however, little of this
potential has been realized.

Bo Ranneby ()
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M.T. Wells and A. SenGupta (eds.), Advances in Directional and Linear Statistics, 201
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Questions related to environmental health, biodiversity, and spatial patterns in
general require new statistical methods. Methods and models must be developed
under so called non-standard conditions — conditions under which many statistical
methods do not work properly but frequently appear in environmental and remote
sensing applications.

In this paper the focus will be on the classification of land use using multispectral
and multitemporal satellite images and some convergence results for nearest neigh-
bour balls (NN-balls). Section 14.2 is devoted to classification methods, including a
brief review of the conventional classification methods, new developments of non-
parametric classification, and the probabilistic classifiers. The reference sample plot
method (today named k-NN) is very popular in natural resource applications. Unfor-
tunately there are no theoretical considerations about the selection of a suitable
metric and the weights. In Sect. 14.3 we will use NN-balls to show how the weights
should be calculated. Some final remarks are given in Sect. 14.4.

14.2 Classification

Supervised classification of remote-sensing images has been widely used as a
powerful means to extract various kinds of information concerning the earth envi-
ronment. The objective of supervised classification in remote sensing is to identify
and partition the pixels comprising the noisy image of an area according to its class,
with the parameters in the model for pixel values estimated from training samples
(ground truths). Usually, the spectral signature is the main aspect of the classes
used to classify the pixels. For multispectral image data acquired by remote sens-
ing devices, one should be aware that they are very complex entities which have
not only spectral attributes (with correlated bands) but also spatial attributes. Proper
utilization of this spatial contextual information, in addition to spectral information,
can improve the classification performance significantly in many applications com-
pared to the conventional noncontextual rules such as discriminant analysis and the
k-nearest neighbour classifier.

A typical problem with the parametric classification methods is the assumption
of (approximative) normality for the feature vector, which is often not fulfilled in
remote sensing applications. For non-normal densities it is nevertheless possible
to use maximum likelihood (ML) classifiers but with other models for the class
distributions. For example, in [18] mixtures of normal distributions are used for
classification of images. Their results indicate a substantial increase in correct clas-
sification rates compared to classification under normal densities. It is worth noting
that mixture models are examples of models when traditional estimation methods
such as ML have a tendency to fail. It has been observed that in remotely sensed
data feature vectors quite frequently possess bimodal or multimodal empirical dis-
tributions. If the training sets are objectively selected empirical distributions of this
kind will become even more common. Therefore general estimation methods, which
give efficient and robust estimates also when traditional methods break down, are
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of fundamental importance for parametric classification. For recent achievements in
this direction we refer to [7, 14, 19].

One way to utilize information from neighbouring pixels is to use the wavelet
transform and denoising techniques. Yu and Ekstrém [23] evaluated various tradi-
tional classifiers by using the wavelet transform on multispectral images, and found
that no matter which type of spatial correlation between pixels the image has and
which kind of conventional classification method one uses, the classification by
using the wavelet transform (specially the wavelet shrinkage) yields a much lower
classification error than that working directly on the original image data, as soon as
we have a class image with some structure in it. It is worth noting that this study is
based on a model for feature vector assuming normality of class distributions.

Concerning the conventional nonparametric classification methods such as the
k-nearest neighbour classifier, its successful combination with the wavelet trans-
form and information theory [15,25] has shown a great potential in remote sensing
applications. The methodology will be described in the following subsections.

We assume that a given pixel from the scene belongs to one of a fixed number of
classes, say, C1, ..., Cg. The proportion of pixels belonging to class C; in the popu-
lation under study is denoted by 7 ;, which is usually unknown. Each pixel gives rise
to certain measurements (e.g., spectral signatures in remote sensing images, or their
various transformations), which form the d-dimensional feature vector X € RY.
Our task is to allocate each pixel into one of the classes in {Cq,...,Ck}, on the
basis of the observed value X = x and/or prior information about the classes.

14.2.1 Conventional Classification Methods

Here follows a short description of three traditional classification methods. See [11,
16] for a comprehensive review of these methods.

14.2.1.1 Parametric Model

In parametric models, the feature vectors from class C; are assumed to be distributed
according to the density p;(x). Then the posterior distribution of the classes after
observing x is p(C;|x) « 1; p(X).

If we assume the probability model in which the observations for class C; are
(d-dimensional) multivariate normal with mean p ;i and covariance matrix X';, the
Bayes rule is to allocate a future observation x to the class that minimises

0; = —2log p(Gj[%) = (x — ;)" 57 (x — ;) + log | Z;| — 2log ;.
This method is known as quadratic discriminant analysis (QDA). When the classes

have a common covariance matrix X', minimizing Q ; is equivalent to maximizing
the linear terms
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Li=2x"Y""'p;, —ul' X7 'pu; +2logm;,

which leads to the linear discriminant analysis (LDA).
In practice, we replace p s Y or ¥, and 7r; with their estimates (e.g., maximum
likelihood) by using a training sample.

14.2.1.2 Nonparametric Model

In nonparametric designs we do not assume any model of the probability distri-
butions. There is a number of nonparametric classifiers based on nonparametric
estimates of the class densities or of the log posterior, such as kernel methods,
orthogonal expansion, projection pursuit, and others. Here we use the simple adap-
tive kernel method which gives a classifier known as the k-nearest neighbour rule.
The k-NN rule is introduced by [6], which is one of the most theoretically elegant
and yet simple classification techniques. It is based on finding the k nearest (usually
in the Euclidean distance) pixels from the training samples (prototypes), and taking
a majority vote amongst the classes of these k samples, or equivalently, estimating
the posterior distributions p(Cj;|x) by the proportions of the classes amongst the k
samples. The nearest neighbour rule (when & is 1) is of special interest in our later
applications.

Nonparametric classification is often associated with the notion “prototype”. A
prototype is a representative point of certain class in the feature space R and is
attached with both field and satellite information. Different classes can have differ-
ent number of prototypes, depending on the property of field data or ground truth
we have. Prototype selection plays a key role in the nonparametric classification.
k-NN is a prototype classifier.

Specifically, assuming the number of prototypes for class C; is n; and n =
Zle nj is the number of all prototypes. For an arbitrary pixel with observed fea-
ture value x, let B(x) denote the hypersphere with centre x and containing exactly
k prototypes and ||B(x)|| the volume of B(x). The number of prototypes in B(x)
from class C; is denoted by k ;. The unconditional probability density of x and the
class-conditional probability density for class C; can be approximated as

k ,
PO e M PN sl (b
respectively. Then the posterior probabilities are obtained as
kj nj
(Gl = L BTEOT 7 _ Ky (14.2)
P) ATB@ k

which implies that the k-NN classification rule assigns the class label independent
of the volume || B(x)||.
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When we are dealing with sparse or rarely occurring objects, however, it is
unrealistic to assume that the number of prototypes for these classes is large so
the approximation above may be violated. Thus there is an obvious risk that such
classes will be underestimated. To overcome this problem alternatives of deriving
the probabilities are needed.

The k-NN classifier uses the whole prototype set of any class to choose the neigh-
borhood, which differs from using the k-nearest neighbour density estimate for each
class. Ties in distances can occur with finite-precision data. One solution to this
problem is to include in the vote all distances equal to the kth largest.

Notice that “nearest” implies a certain metric over the feature space R?. No
matter which metric is used, the k-NN rule is applied in the same way. However,
this metric has to be defined properly because the closeness in the feature space
is no longer so simple as in Euclidean case. This is connected to the similarity in
spatial statistics, which means that the smaller the distance, the higher the similarity
between input data and the prototype.

14.2.2 Nonparametric Classification: New Developments

Very often objects with high environmental values (such as old deciduous forest)
are rare. Remote sensing classification of sparse and rarely occurring objects in
the landscape is complicated and reported classification rates are usually poor. For
several reasons traditional methods will not give satisfactory results:

e The number of field plots in different classes varies too much leading to an
unfavorable treatment of sparsely occurring objects.

e Data editing methods, like the multiedit algorithm [16], often drop whole classes
when applied to moderately sized training sets with more dimensions and classes.

e Noncontextual methods neglect information from neighbouring plots and the
feature vector is not normally distributed.

e Parametric contextual classification methods do utilize information from neigh-
bouring plots but the demands for field data increases, and estimation of some
parameters in the model is unreliable and in many situations impossible.

e The feature vector is not normally distributed and the different class distributions
are often highly overlapping.

To overcome these problems nonparametric approaches have been used. Infor-
mation from neighbouring pixels will be utilized by using the wavelet transform to
denoise the original images by removing small wavelet coefficients and inverting
the wavelet transform. Hereafter a suitable classification algorithm is applied. For
previous work in this direction see [3, 15, 20, 22-26].

The nonparametric classification that we have developed is based on a new
approach for classification of multitemporal satellite data sets, combining multispec-
tral and change detection techniques. The classification method has to be adapted for
handling a mixture of reference data from different sources in order to compensate
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Table 14.1 Definition of forest classes

Proportion deciduous Age
10-39 40-69 70-109 110+
Pine > 70% 1 7 13 19
0-19% Spruce > 70% 2 8 14 20
“Remaining” 3 9 15 21
20-49% 4 10 16 22
50-69% 5 11 17 23
>70% 6 12 18 24

Class 25 = basal area < 1 m*ha™!

for the lack of ground samples from rare classes within the main reference data
source.

As an example, classification of forest land in a study area located in the center
of Sweden is presented. In this application the forest is defined into 24 different
classes with respect to species composition and age, supplemented with the class
“low basal area”, when species composition and age do not have any meaning.
Table 14.1 presents the definition of those 25 forest classes to be used.

The field data from the Swedish National Forest Inventory (NFI) and satellite
images with different dates were used. The dimension of the feature space is 12.

Our classification algorithm is based on the nearest neighbour method. The main
procedures are as follows.

e Define the target function. The probabilities of correct classification for each
class are used, so that each class is of equal importance. Note that it differs from
the overall correct classification, which is often used in various applications.

e Denoise the feature vector. The wavelet shrinkage method based on 2D-wavelet
transform is used to denoise the images. The feature vector consists of compo-
nents that are pixel values from different spectral bands of different temporal
images over both years and seasons.

e Remove outliers from the reference data. When the feature vector has high
dimension and the number of classes is large, new data editing methods are
needed in order to remove outliers due to poor quality of field data, and to find
out the prototypes for each class in the case of nearest neighbour classifiers. This
must be done so that poor quality data is removed at the same time as most of the
natural variations within the different classes are kept. This is achieved by work-
ing with a further subdivision of the classes with respect to other information, as
e.g. basal area, and for the new classes (now 109 classes) suitable techniques for
outlier removal are applied.

e Calculate the information values in the components in the feature vector. Before
the feature vector can be used the components have to be rescaled. The size of
the rescaling factor is a function of the dependence between the studied objects
and the component. Since the classes are defined by more than one variable,
there will not exist any natural order relation between the classes so conven-
tional dependence measures, such as the correlation coefficient, cannot be used.
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Instead measures with the origin in information theory are used. Here we apply
the normed information rate, IRg 5, introduced by [13], which measures the
divergence between the general 2-dimensional distribution and the 2-dimensional
distribution calculated under the assumption about independence and then uses
Shannon’s entropy for the general 2-dimensional distribution to get a measure
between zero (independence) and one (perfect dependence). The definitions are
as follows:

IRe, = 21 (14.3)
where ) )
PE=in=))
PE=i)P(n=))

is the Kullback—Leibler information rate for the class variable & and the feature
component variable 7, and

Iy=> PE=in=j)log

iJ

Hey=—Y PE=in=j)logPE=in=j)
i,j

is the Shannon joint entropy for (&, n).
e Determine a proper metric. We prefer to use the following metric:

d

2
Zwi

i=1

xz(s)q‘xl(t)‘ ) (14.4)

Here x(s) = [x1(s5), x2(8),...,xq(5)], X(®) = [x1(), x2(),...,xq(t)] are two
pixels at location s and ¢, with attribute of the spectral components, d is the
dimension of feature vector, ¢; is the inter-quartile range of the ith feature
component, and the weight

wi = [1 Rclass, feature component (l)]p (14.5)

is determined by the normed information rate of the ith feature component
obtained from (14.3), raised to some power p. And p is determined so the target
function is maximized. Usually p equals to 1.

e Determine prototypes for the classes. Here the information about the occurrence
of different classes in the reference data is used.

¢ Run a nonparametric classification. Here the nearest neighbour classifier is used.

e Declare the quality of classification result by using probability matrices. The
probability matrix is based on the confusion matrix and defined as P = [P;;]
where P;; is the probability that class C; is classified as class C;.

As a result in the example, the probability for correct classification varies
between 30 and 40% for most of the pure classes although it is considerably lower
for some other classes, especially those with different types of mixed forest. The
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Table 14.2 Probabilities of correct classification for C; and Cyy
Class LDA with prior LDA without prior Our method
C 70% 12% 39%
Coy 0% 34% 37%

largest class is C; (younger pine forest) and the smallest is Cy4 (older deciduous
forest). We have compared our classification rates with LDA with or without prior
information. Note that it was impossible to apply QDA here because the field plots
for small classes were too few to be able to estimate the covariances. LDA with prior
information is the most commonly used method. With this method the probability
for correct classification is 0% for 14 out of the 25 classes. The reason is that all
small classes essentially are classified as the larger classes, particularly Cy, which
gets a rate of 70% for correct classification. If we do not use any prior information
then the rates increases for the small classes but drops dramatically (to 12%) for
class C;. A comparison for the largest and smallest classes is given in Table 14.2.

As we want to have high classification accuracy also for small classes they have
to be overestimated (unless the classification is 100% perfect). To overcome this
problem we can calculate matrices for area correction and the precision in the cor-
rection matrices, using Monte Carlo techniques. From this it is possible to obtain
unbiased estimates for the different classes and standard errors of the area estimates.
Resulting estimated areas and standard errors are omitted here.

It is worth to notice that the estimation property is reliable only on the scene
level and it is inappropriate to apply it to the local area or pixel level. To obtain area
estimation of good quality (such as unbiasedness), a new approach is needed, and
we will take it up in the coming subsection.

14.2.3 Probabilistic Classifier

For several applications and a large number of users it is necessary to give pixelwise
information. However, as the distributions for different classes are highly overlap-
ping it is not possible to get satisfactory accuracy at pixel level. It is, therefore,
necessary to introduce a new concept, pixelwise probabilistic classifiers. Instead of
classifying each pixel to a specific class, each pixel is given a probability distribu-
tion describing how likely the different classes are. This new concept has several
important applications: (a) it is possible to give quality declarations at pixel level,
(b) it is possible to derive unbiased area estimates also for small areas and deviating
areas, (c) it is possible to derive cost-efficient monitoring systems.

The concept of pixelwise probabilistic classifiers is closely related to fuzzy clas-
sification; see [2] for different definitions. The drawback with fuzzy classifiers is
that until now the methods are not based on any rigorous theory and the literature is
sparse where the probabilistic classifier definition is used.
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In principle, probabilistic classifiers can be derived both from parametric and
nonparametric classification methods. However, deriving probabilistic classifiers
from parametric methods is a complicated task since for several classes the distri-
butional assumptions are not satisfied. Furthermore, it is of key importance to have
consistent parameter estimates. To avoid the problem of misspecification, nonpara-
metric approaches are preferred. The probabilistic classifier derived here is based on
the k-NN method with k = 1, defined in Sect. 14.2.1.2.

Articles on fuzzy classification and k-NN are available, see e.g. [1, 10], but they
have a different scope. To be useful for area estimation and quality assessment at
pixel level the probabilities must be extracted from proper probability distributions
for the different classes. When the classifier uses distances in feature space to derive
the probabilities it is intuitively obvious that the probabilities will be inversely pro-
portional to the distances raised to some power. However, there is only one value
that is correct and that value depends on the dimension of the feature vector. Actu-
ally, by using the nonparametric density estimation [12], the density for class C; atx
will be inversely proportional to the volume of the nearest neighbour ball with cen-
ter x. Note that the volume is proportional to the radius, based on the metric defined
in (14.4), raised to the dimension of the feature space. The derivation is similar as
for (14.1) and (14.2), but now using the NN-balls in the feature space.

As a theoretical background, we introduce a proposition based on [14] before the
derivation. Let B(x,r) = {y : |[x —y| < r} denote the ball of radius r with center

at x in the feature space. Then the volume of the ball B(x,r) is || B(x,r)| = cqr?
where cqg = 7%/2/I'(d /2 + 1).
Let X1,X>,..., X, be a sequence of i.i.d. d-dimensional random vectors with

the common density function g(x) and define the nearest neighbour distance to the
point X; by
Ry (i) = min [X; — X;|.
J#i

Then B(X;, R,(i)) is the so-called NN-ball of X;.

Proposition 14.1. Let X1,X5,...,X,, be i.i.d. random vectors with the common
density function g(x) and

Y (i) = n ||B(X;, Ru(i))|| = nca R4 (i).

Then b
X;, Y,(0) — (X,Y), asn— oo, (14.6)

where X has density g(X) and Y given X = X has an exponential distribution with
parameter g(X).

Note that (X;,Y,(i)),i = 1,2,...,n are exchangeable and asymptotically
independent, and the joint probability density of (X, Y) is g2(x) exp{—yg(x)}.
Furthermore,

E[Y,()X; =x] > E[Y[X =x] = 1/g(x).
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Let now

R, (x, C;) = the minimum distance in the feature space (using the metric (14.4))
from x to the prototypes of class C;. 14.7)

The conditional probability density for class C; can be estimated by

1
pj(x) = . (14.8)
! nj | Bx. Ru(x.C)))|
Then the posterior probabilities are obtained as
R m;pj(x)
PO =
Zi:l i pi (X)
T Z T
n; HB(X, R,,(X,C-))” Pl | B(x, R, (x, C;))|
w; _n]/n Z
- nmxmuC»n _JMxm@cm
= n " (X’ CJ) .
Yini Ry (x.Ch)
For each pixel with location s and feature vector x(s) = [x1(s), x2(s), ..., xg ()],

we assign the following proper probability to class C;

: R4 (x(s), C))
I(s) = L . 14.9
re Y RyA(x(s), Cr) (142

Thus, the probability vector at pixel s is obtained as p(s) = [p'(s). p?(s).....
K
PG

14.2.4 Applications of the Probabilistic Classifier

The probabilistic classifiers can be used in different ways. For instance, it can be
used to measure uncertainty at pixel level, or to obtain reliable area estimates locally.
It can also be applied to correct the erroneous mask of maps (see comments in the
last section).

e Quality evaluation of traditional classification
A traditional classification is obtained by choosing the class with the highest
probability for each pixel. With the probabilistic classifier, it is possible to give
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quality declaration at pixel level. The pixelwise vectors of probabilities (14.9) can
be used to judge how reliable a traditional classification is and to derive measures
of the uncertainty (entropy) for the individual pixels. Based on the probability
vector, the entropy at pixel s is defined as

K
- Z P’ (s) log p’ (s). (14.10)
j=1

e Monitoring systems
In application, in addition to the classified map a raster map expressing the qual-
ity (entropy defined in (14.10)) for each pixel is produced. Areas where the
precision is unsatisfactory will be identified. In these areas it is possible to take
additional field plots and perform an improved classification. Here it is important
that the classification method can handle different types of reference data. The
final product will be two maps, one showing the pixel wise classification and
the other giving the classification accuracy at pixel level. Figure 14.1 illustrates
our concept of probabilistic classifiers operated on a small site of size 16 x 16
pixels. The area consists of mixed forest, which implies difficulty to classify.

E:

xample of entropy and probability calculation

%
81 82 83 84 85 8 87 8 89 90 91 92 93 94 95 96
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egend - entropy
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[ 0.75-1
1-1.25
1.25-1.5
1.56-1.75
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. E2-225
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w1 Field
el o ]
< investigations in !

i selected areas L

Fig. 14.1 The concept of probabilistic classifiers. Test area with mixed forest, eight classes. Field
plots can be added in areas where entropy is high (i.e., quality at pixel level is unsatisfactory) to
produce additional reference data as input to a renewed (improved) classification
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Darkest colored pixels indicate higher entropy values or unsatisfactory quality.
The diagram on top right shows the probability distributions of 16 pixels (one
row) picked from the entropy map on the left side. For a given pixel, the more
even distributed probabilities among classes, the high entropy. If one class domi-
nates the probability vector (i.e., having high probability), then the pixel has very
low entropy and is reliably classified.

Area estimation

Although it is possible to obtain satisfactory area estimates on scene level using
area correction matrices derived from the confusion matrices as we discussed in
Sect. 14.2.2, the precision is not good enough for smaller areas or on pixel level.
The pixelwise vectors of probabilities can be used to get unbiased area estimates
also for quite small areas and for deviating areas, such as potential areas for
nature reserves. The area estimates is obtained simply by summation of the “class
probabilities” over the selected subregion. It is extremely important that proper
probability distributions allowing frequency interpretation are derived; otherwise
misleading results are obtained.

Concerning the quality assessment related to our new area estimates, resam-
pling and subsampling methods will be considered. The advantage of such
methods is that statistical inference can be done without knowledge of the under-
lying spatial dependence mechanism and marginal distributions that generated
the data. Furthermore, for the user no explicit theoretical derivation is necessary
but instead intensive computing. Most of the proposed methods in the literature
assume stationary data. However, when modeling real-life data, the hypothesis
of stationarity often must be rejected. Especially for nonstationary spatial data,
there are very few available methods to do inference. Sherman [17] has provided
a short discussion on subsampling of spatial lattice data in the context of regres-
sion models with stationary errors, and later [4, 5] has proposed resampling and
subsampling methods for nonstationary spatial lattice data. The nonparametric
variance estimator proposed by them can be used to estimate not only the vari-
ance of the area estimates derived from the probabilistic classification, but also
the variance of estimators for continuous variables (such as wood volume) with
respect to (estimated) area of interest (e.g., forest).

14.3 k-NN Estimation in Natural Resources

Starting around 1990 with articles by [9, 21], k-NN techniques have gained pop-
ularity for natural resources applications. Kilkki and Pivinen [9] used the term
“reference sample plot method”. Later the term “k-NN method” was established
although the method differs from the k-NN method introduced by [6]. Katila and
Tomppo [8] provided a survey of the early k-NN (reference sample plot method)
literature for estimating forest attributes. For prediction we have observations of
the response variable Y for a subset of pixels and observations of covariates X
(d-dimensional feature vector) for all pixels. The k-NN prediction for the i th pixel is
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-1
k k
Z Wij Z WijYij»
j=1 j=1

where {y;;, j = 1,2,...,k} is the set of observations closest to the ith pixel with
respect to a metric in the feature space, and w;; is the weight assigned to the jth
nearest neighbour observation. Common selections for the metric are Mahalanobis
distance or Euclidean distance. Usually the weights are chosen to be inversely pro-
portional to the distance raised to the power of 1 or 2, regardless of the dimension
of the feature space.

Theoretical considerations about the selection of the weights and a suitable met-
ric are lacking. For selection of a suitable metric some of the ideas described in
Sect. 14.2.2 will be useful. In this section we will use NN-balls to show how the
weights should be calculated.

For the reference set (e.g., satellite data) we have information both for the
response variable Y and the feature vector X. For a pixel with value x, the best pre-
dictor of Y, given squared loss function, is the conditional expectation E[Y |X = x].
It can be written as

E[Y|X =] = [ (v Ix)dy = / y%w (14.11)

The support of Y is partitioned into sets C;, j = 1,..., K. Then

Ko
EYX=x]=% — d
[Y[X =x] ;p(x)/ciyp(xw)p(y)y
K
1 yp(y)
=N ___pyec) | 22 dy.
Lo 0w e e

If the partition {{_J C;} is fine enough then p(x|y) is almost constant for all y € C;,
and denoted by p(x|C;). This gives us

K

1
B == 3Pt € Gy [ s
J=1 i j
= Z (X)P<Y € C)pXIC)1;. (14.12)

Here 1 ; is the expected value of the response variable for class C; and p(x|C;) is
the density of the covariates for class C;.

Suppose now that for the class C; we have n; reference points and let n =
Zle nj, the total number of reference points. Suppose that a suitable metric in
the feature space, such as (14.4), has been chosen. With respect to that metric the
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distance from the pixel with covariate value x to the nearest pixel in the reference
set belonging to class C; is denoted, as in (14.7), by R, (x, C;). As we have seen
before the volume of the nearest ball converges in distribution to an exponential
distribution,

nj; ”B(X, Rn(X, Cj)) ” = I’ledRZ(X, Cj) 2) Zj,

where Z; is exponential distributed with mean 1/ p(x|C;). Furthermore,

1

EnjcqRI(x.C))] — E[Z;] = PR
]

To obtain a predictor y(x) of the pixel with covariate value x we substitute each
component in the best predictor E[Y |X = x| (14.12) with a reasonable estimate.

Thus,
K

. 1
yx) = Z S0 ¥ € CAHICHA,. (14.13)
If the reference set is chosen properly, then
nj
— — P(Y € Cj).
n
The mean pt; has to be estimated by the observed value y ; Vi and the density p(x|C; )

will be estimated by [1 ;¢4 Rd (x,C;)]™1, and p(x) by Z [njcdR (x,Cp)]!
Thus

_ln

yx) = p(x) Zyj—[n]cdR x G

K
=Y Rx.C) Zy, R;4(x.C)). (14.14)

Consequently, the weights in k-NN estimation shall be inversely proportional to the
distance raised to the power of d, the dimension of the covariate (feature) space,
and not to 1 or 2 which are standard choices in the k-NN (reference sample plot
method) literature.

14.4 Final Remarks

Remote sensing classification of sparse and rarely occurring objects in the land-
scape is complicated. An approach, based on nonparametric statistical methods,
has been developed to overcome the problems arisen with traditional classification
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methods. It utilises information from multitemporal and multispectral satellite data
and change detection techniques. The classification method is also adapted for han-
dling a mixture of reference data from different sources in order to compensate for
the lack of ground samples from rare classes within the main reference data source.
The new algorithm is derived in order to optimize the average probability for correct
classification, i.e., each class is equally important.

As we have seen the probabilistic classifiers are useful for measuring uncertainty
at pixel level and obtaining reliable area estimates locally. It can also be applied
to correct the masks of maps. The masks from the topographic maps are usually
used to define different land use of vegetation, e.g., forest land, peat land, or agri-
cultural land. Then the classification is performed within the mask. It has, however,
been found that the errors in masks can be significant, and thus development of bet-
ter masks is needed. Otherwise we know a priori that the classification accuracy
will not be satisfactory. An application of the probabilistic classifiers to obtain new
masks for forest land and peatland has been conducted successfully in other studies
of the authors. The idea is as follows: (a) use the probabilistic classifier together
with information from digital elevation model (wetness index) and neighbouring
pixels to get the probability vector for classes (both forest and peatland) at pixel
level and consider it as a prior probability distribution for each pixel; (b) apply the
probabilistic classifier by using remotely sensed data to get the probability vector
for classes at pixel level; (c) join (a) with (b) to get the a posterior probability vector
for each pixel; (d) aggregate the classes to two categories: Forest and Peatland; (e)
calculate entropies for each pixel and determine the threshold value according to
the entropies so that pixels belong to one of the following zones: Forest, Peatland,
or Transition Zone; (f) recalculate the probability vectors for pixels in Forest and
Peatland zones. In such a way, two masks with higher quality are obtained for forest
and peatland, respectively. A third one, the Transition Zone, is given for pixels with
a certain degree of uncertainty.
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Chapter 15
Probabilistic Recurrence Relations

M. Bhaskara Rao, S. Kasala, and H. Zhang

Abstract A sampling of discrete probability problems, some of them coming from
consulting work, is presented. We demonstrate how a probabilistic recurrence rela-
tion arises from the pit of the problem and present ways and means of solving the
recurrence relation.

15.1 Introduction

Recurrence relations crop up frequently in mathematical research. The basic set-
up is as follows. A sequence X,,n > 0 of numbers is said to satisfy a recurrence
relation of order k > 1 if

Xp=a1Xna+aXp o+ +arXnk +bp.n>k

for some fixed constants aq,as,--- ,ag, and sequence b,,n > k. The goal is to
find an explicit mathematical expression for the nth term X,. In order to find an
expression, we need the values of Xo, X1, , Xr—1. These values are collectively
called ‘boundary conditions’. The mathematical form, of course, depends on the
‘boundary conditions’. If it is not possible to find an explicit mathematical expres-
sion for the nth term of the sequence, one could investigate the asymptotic behavior
of the sequence. If the sequence b,,n > k is a constant, i.e., b, = b foralln > k,
the recurrence relation is said to be homogeneous. If the relation is homogeneous,
one could use ‘difference equations’ methodology to find an explicit expression for
the nth term of the sequence. If the relation is not homogeneous, one has to devise
some ‘ad hoc’ method to solve the recurrence relation.
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A classical example of a recurrence relation is the one satisfied by the Fibonacci
sequence X,,n > 0,ie., X, = X,—1 + Xn—2,n > 2 with boundary conditions
Xo = 0and X; = 1. This is a homogeneous recurrence relation of order 2. The nth
term has an explicit expression:

_ 9" -1 —-9)"

X, = n >0,

N

where ¢ = @ is the so-called Golden Ratio.

In this article, we focus on recurrence relations that arise from probabilistic
investigations. Probabilistic recurrence relations are of two kinds. The sequence
Xn,n > 0 could be a sequence of probabilities satisfying a recurrence relation
of order k for some k > 1. Solving such recurrence relations falls into the realm
of mathematics. On the other hand, the sequence X,,n > 0 could be a sequence of
random variables satisfying a recurrence relation of the type

d
Xn = f(Xpn—1, Xn—2,.... Xpn—r), n=>k,

where the symbol 2 tmeans ‘equal in distribution’ and the distribution of X, is the
same as the distribution of f(X,—1, Xn—2,..., Xy—). The function f(-) is fixed
and known. Typically, the function f(-) is of the form

f(Xn—h Xn2,..., Xn—k) =a1Xp-1+ax Xy o+ -+ apr Xy + Yy,

where ay,as,...,ay are constants and Y, is a sequence of random variables inde-
pendent of the sequence X,,n > 0. The goal is to determine the distribution and
properties of X,,. In order to determine the distribution and properties of X, such as
mean and variance, we ought to have a knowledge of the distribution of the initial
random variables Xg, X1, ..., Xx_1, and also that of Y;,,n > k.

The probabilistic recurrence relation outlined above looks like an Autoregressive
model encountered in Time Series Analysis but the random variables involved have
special distributions. In our ‘modus operandi’ the random variables have discrete
distributions. Further, we do discuss situations in which the recurrence relations are
not exactly the type outlined above but in spirit they are similar.

In this article, we present a number of examples from discrete probability which
are unified by the presence of a probabilistic recurrence relation. Some of these
problems came from our consulting work. These are listed below:

Dermal Patch Problem
Patterns in Coin Tossing
Chemical Bonding Problem
Yell Game

Noodles Problem

Nk e =
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These examples can be used in a class-room setting to demonstrate how proba-
bilistic recurrence relations arise naturally in some probabilistic investigations and
exhort students to find ways to solve such recurrence relations.

15.2 An Illustrative Example

Let X1, X5, -+ be a sequence of independent identically distributed Bernoulli ran-
dom variables with Pr(X; = 1) = p =1—Pr(X; = 0) with0 < p < 1 fixed.
What is the distribution of S, = X; + X5 + --- + X,,? There are many ways this
problem can be solved. We can solve this problem by building a probabilistic recur-
rence relation. Note that S, = S,—; + X,. We postulate the induction hypothesis
that S, ~ Binomial(n, p) foralln > 1,i.e., Pr(S, =r) = ('r')p’(l -p)"r=
0,1,---,n. The result is true for n = 1. Assume that the result is true for all k < n.
We will show that the result is true also for k = n + 1. Observe that S, 4+, = 0 if
and only if S, = 0 and X,,+; = 0. In view of independence,

Pr(Sys1=0)=Pr(S, =0)-Pr(X,41 =0) = (1 - p)"t1

Let 1 <r < n.Observe that S, = r if and only if S,, = r and X,,4+; = O or
Sp =r —1land X,,4+1 = 1. In view of independence,

Pr(Sy+1=r)=Pr(S, =r)-Pr(Xy+1 =0 +Pr(S, =r—1)-Pr(X,+1 =1)

B (lj)”r(l —-p)" - (=p)+ (, " 1)19"‘(1 —p) T
()L o
r r—1
1
) <+ )”r“—pr’—rﬂ

Finally, S,,+1 = n + 1 if and only if S,, = n and X,,+; = 1. Consequently,
Pr(Sps1=n+1)=Pr(Sy =n)-Pr(Xps1 =1)=p"-p=p"tL

Thus S, +1 ~ Binomial(n + 1, p).

15.3 Dermal Patch Problem

A dermal patch has been created to release doses incrementally over a period of
10days into an organism to which the patch has been attached. The goal of the
doses is to reach ten receptors, currently dormant, inside the organism, and make
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them active. On Day 1, the patch releases one dose, which latches onto one receptor
and makes it active. On Day 2, the patch releases two doses, which latch onto two
receptors, one dose per receptor. If the receptor is already active, the new dose makes
it inactive. If the receptor is not active, the new dose makes it active. On Day 3, the
patch releases three doses of medicine, which latch onto three receptors, one dose
per receptor. The active or inactive status of the receptor which received a dose
follows the same paradigm outlined above. This will continue until the tenth day,
on which ten doses are released, which latch onto all the ten receptors, one dose per
receptor. The question, posed by a pharmaceutical company, is to determine how
many receptors are expected to be active at the end of the tenth day.

We have generalized and synthesized the problem. We have christened the
generalization as the ‘Light Bulb’ problem.

In the light bulb problem, we assume that there are n > 1 light bulbs, serially
numbered from 1 to n, each one attached to a toggle switch. If the bulb is off and
its switch is pressed, the bulb lights up. If the bulb is on and its switch is pressed,
then the bulb is off. Initially all bulbs are set off. On Day i(= 1,2,...,n) exactly
i of the n switches are randomly pressed. The question raised is to determine the
expected number of light bulbs on at the end of the nth day. The problem was first
considered in [8].

Let X, be the number of light bulbs on at the end of day n. Write

Xn=Xum + Xuz + -+ Xy,

where X,; = 1,if the ith bulb is on at the end of Day n, and = 0 otherwise. Note that
Xn1, Xn2,- -+, Xnn are exchangeable. It suffices to calculate pi") =Pr(X,; =1).
This would give E(X,) = np™. Let

pir) = Probability that the bulb attached to Switch 1 is on at the end of Day r.
Then the light bulb attached to Switch 1 lights up at the end of Day r if and only if
(1) the light bulb was on at the end of Day (r — 1) and Switch 1 was not pressed on
the rth day; or (2) the light bulb was off at the end of Day (» — 1) and Switch 1 was

not pressed on the rth day. We then come up with the following recurrence relation,
a non-homogeneous difference equation of order 1,

r _ r _ r 2r _
p = (=) S l))=—+(1——)pir Vor=23 .,
n n n n

r)_1

with boundary condition pgl) = 1/n. To solve this equation, letting qﬁ” =p =3

r =1,2,---,n, we have the following recurrence relation

0 2r\ -1
=(1-= i
q1 ( n )‘11
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forr = 2,3,.--, n with boundary condition qgl) = % — % Thus, we obtain
o 1 (1 1\{ 2i 1 d 2i
W=+ (30 7) :z(l‘n(l‘z))’
i=2 i=1
forr =1,2,---,n. Therefore,

E(Xn)=%(1—ﬁ(1—%)).

i=1

Note that £X,, = g if n is even, and ~ % if n is odd. The idea of recurrence relation
has been extensively exploited in [8] to calculate the variance, covariance and the
distribution of Xj,.

15.4 Patterns in Coin Tossing

If we toss a fair coin four times, what are the chances of getting the pattern HHHH?
HHHT? They are the same. Let us pose the problem differently. Keep tossing the
coin until we get the pattern HHHH. How many tosses are needed to get the pat-
tern HHHH? How many tosses are needed to get the pattern HHHT? It is harder to
get the pattern HHHH than the pattern HHHT in the sense that the expected num-
ber of tosses required to get the pattern HHHH is greater than the corresponding
expectation for HHHT.

Questions on expected number of tosses needed for any pattern of outcomes to
appear for the first time have been solved elegantly using martingale arguments
in [6] and [5]. Their work was further extended in [7]. In this section, we focus
on the distribution of the number of tosses required. We show how probabilistic
recurrence relations help solve the problem and provide a rich source and variety of
number sequences.

Let Ty be the number of tosses required to get heads for the first time. It is an
elementary exercise that the distribution of Ty is geometric, i.e.,

Tu: 1 2 3
Probability: 12 (1/2)2  (1/2)3

with E(Ty) = 2.
Let Ty be the number of tosses required to get the pattern HH. It is not hard to
show that its distribution is given by

Probability: (1/2)2 (1/2)3 2(1/2)* 3(1/2)° 5(1/2)°
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with E(Tyg) = 6. A surprising feature about the distribution is that the coeffi-
cients of power of 1/2 form the Fibonacci sequence! The distribution of Ty can be
derived from that of Ty. The key idea is a probabilistic recurrence relation.

Suppose that we are observing Ty. Look at the (T + 1)® toss. It could be either
H with probability 1/2 or T with probability 1/2. If it is H, we have the pattern
HH occurring for the first time and Tyg = 1 + Ty. If it is T, we need to start
from scratch beyond the (Ty + 1)* toss to look for the pattern HH. Consequently,
Tun = 1+ Tu+Tijy. where Ty is an independent copy of Tuy. These deliberations
lead to the following probabilistic recurrence relation

1+ Ty with probability 1/2,

Toy =
i { 1 + Ty + Ty with probability 1/2.

It is easy to exploit this recurrence relation to get E Tyy.
1 . 1
ETgg = (1 4+ ETy) - 3 + (1 + ETyn + ETyy) - 3
=3+ ! ET;
= ) HH,

implying that ETyy = 6. This relation can be exploited to calculate variance of Tyy
and distribution of Tyy. As for the distribution of Ty, note that

Tyg =2 ifandonlyif Ty =1.

Consequently,
P(Th 2) ! Pr(T; 1) hy’
= = — r = = — .
HH 3 H >
Also,
Tyn =3 ifandonlyif Ty = 2.
Hence,
P(Tyg =3) = ! Pr(Ty=2) = 1y’
HH = 2) = {5 rn = 2) = 7] -
Now
Tyu =4 ifandonlyif Ty =3 or
THH =1 and TI:H =12.
Therefore,

-(3) () 6) () =2()
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One can proceed inductively to lay out the entire distribution of Tyy. It should be
pointed out that there are different ways to develop a recurrence relation for Tyy.
Following the same trail, we have obtained the distribution of Tyyy.

Tyun: 3 4 5 6 7 8 9
Probability: (1/2)® (1/2)* 2(1/2)5 4(1/2)5 7(1/2)7 13(1/2)8 24(1/2)° ---

A serendipitous discovery is that the coefficients of powers of ( %) in the distri-
bution constitute a tribonacci sequence, i.e.,

fi=1Lfa=1 =2 fa=4 fs=7, fe =13,
f7 = 24,"' ,fn = fn—l +fn—2+fn—37

etc. In general, one can show that the coefficients of powers of (%) in the distribution
of Ty ... g constitute a n-bonacci sequence. Further,
——

E(Tgy...g)=2"""-2.
N’

Using similar ideas, we have obtained the distribution of Tyr.

Tyr: 2 3 4 5
Probability:  (1/2)2  2(1/2)*  3(1/2)*  4(1/2)5

with E(Tyr) = 4. Note that the coefficients of powers of (%) in the distribution
constitute the entire sequence of natural numbers.

A very rich family of distributions on {0, 1,2, 3,--- , } can be generated by look-
ing at the distributions of Tpaem for a variety of patterns. Further, by looking at
the coefficients of powers of (%) in the distributions, we will have a rich variety
of positive integer sequences. In [4], expectations for patterns of length 3 are given
using Markov Chain ideas. Here is a partial list of expectations.

Pattern Expectation

H 2
HH 6
HT 4

HHH 14
HHT 8

HTH 10
THH 8

One can provide a general formula for E(Tpauen) for any given ‘Pattern’ of heads
and tails. If the ‘Pattern’ is of length n, then
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E(Tpattem) = 2n + bl ° 2n—1 + bZ * 2n—2 + b + bn—l ° 2’

for some by, by, -+, by—1 in {0, 1}. The following algorithm gives the coefficients.

Letaja; - --ay be a pattern of length n.

Stage 0:  Write the pattern as follows.

ayas---dy

aias---dp

There is a perfect alignment.

Stage 1: Slide the second pattern one position to the right.
aijdads---dpy
apdaz---dp—14dn

If there is a perfect match in the common positions of both the patterns, b; = 1,

otherwise, b; = 0.
Stage 2:  Slide the second pattern two positions to the right.

aijdadsdyg - -dy
apdz - --dp—2dn—1dn

If there is a perfect match in the common positions of both the patterns, b, = 1,

otherwise, b, = 0, andsoon .. ..
As an illustration, look at the pattern HHTTHH.

Stage 0:  Write the pattern as follows.

HHTTHH
HHTTHH

There is a perfect alignment.
Stage 1: Slide the second pattern one position to the right.

H HTTHH
HHTTHH
b1 =0.
Stage 2:  Slide the pattern two positions to the right.
HH TTHH
HHTTHH

by

Il
o
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Stage 3: Slide the pattern three positions to the right.

HHT THH
HHTTHH
b3 =0
Stage 4:  Slide the pattern four positions to the right.
HHTT HH
HHTTHH
by =1
Stage 5:  Slide the pattern five positions to the right.
HHTTH H
HHTTHH
bs =1
Consequently,

E(Tunrran) = 2% + 2% +2' = 70.

15.5 Chemical Bonding Problem

A certain system has ten molecules in some hierarchical order from 1 to 10, say. A
catalyst is added to the system and the resultant reaction lines up the molecules. One
can read the hierarchies of the molecules from left to right. If there are molecules in
the line-up in consecutive increasing hierarchical order, then these molecules merge
to become one molecule. A new hierarchical order sets in among the molecules that
are left over including the merged ones. Again, a catalyst is added to the system and
the whole process of merging renews again. The process continues until we get a
single molecule. The question of interest is how many times catalysts are expected
to be added until we get a single molecule.

We have generalized and synthesized the problem. We relabel the problem as
‘Card Shuffling Problem’. (A related problem has been presented in [2]).

In the card shuffling problem, we assume that n cards, serially numbered from
1 to n, are shuffled thoroughly and spread out in a line on a table facing up. The
cards were then read from left to right, and those cards with consecutive numbers
in increasing order are merged. After the mergers, the cards are renumbered serially
from 1 onwards to whatever number of cards available now. Cards are shuffled thor-
oughly again and laid out on a table in a line facing up, etc. The process continues
until only one card is left. See [9] for more details.
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Let X, be the number of shuffles needed so that there is only one card left at
the end. The principal questions are: E(X})? Var(X,)? Distribution of X,,? We will
start with a special case n = 2 to illustrate our approach to the problem, and then
extend the idea to the general case.

Assume n = 2, let )22 be an independent copy of X5. X, takes values {1, 2,
3, ---}. If the first shuffle ends up with the permutation 1 2 (with probability 1/2),
only one shuffle is needed. However, if the first shuffle ends with the permutation 2 1
(with probability 1/2), then the cards are needed to be shuffled again. The number
of shuffles needed to have one card left is X,. In summary, we have the following.

Possible permutations: ~ Probability =~ Value of X,
{12} 1/2 1
21 1/2 14 X,

In other words,

X, — 1 with prob. 1/2, corresponding to the shuffle result {1 2},
2711+ X, with prob. 1/2, corresponding to the shuffle result {2 1},

This is a probabilistic recurrence relation. It can be exploited to calculate E(X>).
For example

E(G) = 16 (1/2) 4 B+ K) % (1/2) = 1+ SE(Xa),

implying that E(X,) = 2.

Before moving to the general case, we follow the notation from [2, p. 168], by
letting T, s be the number of permutations of {1,2,---,n} such that in each per-
mutation there are s pairs of consecutive increasing integers in the permutation. For
n < 8, Ty is given by Table 1 on the same page. We further let A(n, k), k =
1,2,---, n be the number of permutations of {1,2,---,n} in which k cards are left
after merging from n cards. It is shown in [9] that A(n,k) = T(n,n — k) and
Y ko1 A(n k) = n!.

Suppose that there are n cards initially. Let X be an independent copy of X,
k = 2,3,---,n. If the first shuffle ends up with the permutation 12 --- n (with
probability A(n, 1)/n! = 1/n!), only one shuffle is needed. In general, if there are
k cards left (with probability A(n,k)/n!, k = 2,3, ---) after the first shuffle and all
possible mergers, then the k cards are needed to be shuffled again and the number
of shuffles needed to have one card left is X k- Therefore, we have the following
recurrence relation.

1 with probability A(n, 1)/n!,
Y. — 1 + X, with probability A(n,2)/n!
n =

1 + X, with probability A(n,n)/n!
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Hence, we have

A(:;Z)E(Xz) P An,n — I)E(Xn—l) L A(n,n)

E(X,) =1+ E(Xn).

n! n!

and so

n—1
E(X) = (1 £y A(Z;k)E(Xk)) /(1-42).
k=2 ' '

The following table gives E(X,) when n < 10. More results can be found in [9].

n 2 3 4 5 6 7 8 9 10
E(X,) 2 333 461 584 7.02 817 930 1042 1140

15.6 Yell Game

This game is played by many improved comedy troupes for encouraging coopera-
tion between performers. See [1] for details. The game is played by N individuals
who stand in a circle, initially looking down at the floor. On a signal, all play-
ers simultaneously lift their gaze from the floor and each player fixes his/her gaze
exactly at one other player in the circle. If any two players make eye contact with
each other, they yell at each other and they are out of the game. The game continues
with players who remain. In [1], the focus was on calculating probability of silence.
In this section, we focus on determining the distribution of the number of people
left over at each turn of the game.

Let n>2. Let XO(") =n = the number of players at the start of the game. Let
X IE") = the number of players at the end of kth turn of the game. If n is even,
the possible values of X,E") are 0,2,---,n. If n is odd, the possible values of
X,En) are 1,3,.--,n. Let us look at some special cases. Suppose n =2. Then

Xéz) =2, Xl(z) =0, X2(2) =0,--- etc. Suppose n = 3. The distribution of XI(B) is
given by

x®: 1 3
Probability 3/4 1/4

It is clear that X 153), k > 0 constitutes a Markov Chain with state space {1, 3}
and transition probability matrix

F= (3}4 1(/)4)'
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Using Markov Chain ideas or a recurrence relation approach, one can show that the
distribution of X 153) is

@),
xP: 1 3
1

Probability - L

I

with E(X ;3)) =1+ %k. Let us look at the case n = 4. The distribution of X 1(4) is
given by

x®. 0 2 4
Probability 1/27 8/27 18/27

and E(X 1(4)) = 88/27. The sequence X 154), k > 0 constitutes a Markov Chain with
state space {0, 2, 4} and transition probability matrix

1 0 0
P=| 1 0o o0
1/27 8/27 18/27

Using Markov Chain ideas or a recurrence relation approach, one can show that the
distribution of X ]§4) is

x®: 0 2 4
. k. k. k.
Probability 11— g T JS’ 32k +42 32k +92
with E(X 154)) = 2];:12” . We do not have a general formula for the distribution of

X IE") for any n. One interesting problem is to find an expression for the probability
of ‘silence’ for any 7. More precisely, let

pn =Pr(X l(n) = n) = Pr(all players stay in the game at the end of the first turn)
= Pr(No yells).

It will be interesting to obtain an asymptotic formula for p,,.

15.7 Noodles Problem

15.7.1 Introduction

There are n strings. Each string has two ends. Out of 2n ends two ends are selected
at random and tied. If the two ends come from the same string, we will have a loop
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right away. Otherwise, we will have (n—1) strings with 2(n—1) ends. We now select
two ends at random from these 2(n — 1) strings. The selection will continue until
the last two ends available are tied. Let X,, be the total number of loops that will
form. The possible values of X, are 1,2,---,n. The objective of this section is to
determine the distribution of X, its expected value, the variance, and the asymptotic
normality.

15.7.2 Expected Value

The random variables X, and X, have a connection. If the initial selection of
two ends come from the same string, we will have a loop right away and X, =
1 + X,,—1. On the other hand, if the initial selection of two ends come from two
different strings, then X, = X,_;. The probability that two ends come from the
same string is given by 1/(2n — 1). The complementary event has the probability of
2(n — 1)/(2n — 1). Formally, the relationship between the random variables can be
written as

d {14+ X,—1 withprob. 1/(2n — 1),
Xn = 15.1
" Xn—1 with prob. 2(n — 1)/(2n — 1). ( )

This fundamental recurrence relation in distribution can provide formulas for
E(Xy) and Var(Xy). In addition, the distribution of X, is derivable. First, let us
work with the expected value. Observe that

1 2(n—1
E(Xn) = Ed+ X+ 2=V ex, )
2n —1 2n —1
1
= E(X,— =2.,3,---.
2n—1+ ( n 1)»” , D,

The boundary conditionis £(X;) = 1. The formula for the expected value can be
set out directly from the recurrence relation between two consecutive expectations.

E(X)—1+1+1+ + !
Y305 2n—1
Note that the expected value diverges to oo, rather slowly, at the pace of Inn. An
approximation for the expected value is given by

Inn 1
where y ~ 0.5772 is the Euler’s constant. This approximation is good with agree-
ment between the exact value and the approximation taking place in the first two
decimal places for n equal to as low as 15.
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15.7.3 Variance

The recurrence relation between the random variables X, and X,,—; can be exploited
to get a recurrence relation between Var(X,) and Var(X,—1). However, we present
another relation between X, and X,—; by introducing a Bernoulli random variable.
In the context of n strings, let

7 = 1 if the randomly chosen ends come from the same string, (15.2)
"7 | 0 otherwise. '

The probability distribution of I, is given by

P(I, =1)=

=1-P(I, =0).
2n—1 (I =0)

The distributional recurrence relation (15.1) is equivalent to

Xo L Xoot + 1 (15.3)

with X, _; and [, independently distributed. Consequently, in view of independence
of X,,_1 and [,

2(n—1
Var(X,) = Var(X,—1) + Var(ly) = Var(X,—1) + —(2(n 1))2,11 =2,3,---
n —

The boundary condition is given by Var(X;) = 0. The formula for the variance
works out to be

Var(X,) lhipli 1 LI I
ar e - - j— J— J— S .
g 35 2n—1 32 1 52 (2n —1)2

The variance also diverges to oo, rather slowly, at the pace of In . An approximation

to the variance is given by

Var(X,) = 2 {1n2 4 ) 3
ar =—+1In —y -
n = 2" 46

15.7.4 Distribution

The recurrence relation (15.1) and (15.3) can be used for obtaining the distribution
of X,, from that of X,,_;. Let the distribution of X,,—; be given by
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)(n—l 1 2 cee n __1
Probability Pn—1,1 Pn—1,2 e Pn—1,n—1-

Let us denote the probability distribution of X,—; by the (n — 1)-tuple: (pn—1,1,
Pn—12.""* s Pn—1,n—1). From (15.2), following the same notation, the probability
distribution of X, is given by

(pn,h Pn2,:°°, pn,n) = m—1 * (0, Pn—-1,1,Pn—1,2,""", pn—l,n—l)
2n —2

+2n—1

* (Pn—1,1, Pn—=1,2,""* » Pn—1,n—1,0).

The distribution of X, is a convex combination of two probability n-tuples created
from the distribution of X, _1. From this, it follows that, forn = 2,3, ---,

2n —2 2n—-2)2n—4)---(H(2)
Pn1 =5 Pr11 = 2n—1)2n—=3)---(503)’
1 2n—2 .
Pn,j = mpn—l,j—1 + mp”_l’j’] =2,3,---,n—1;
_ 1 _ 1
Pnn = o — 1pn—l,n—l = 2n —1)(2n _3)“.(5)(3).

The distribution of X, for selected values of n is tabulated below.

The distribution of X},
n 1 2 3 4 5 6
1 1 0 0 0 0 0
2 2 3 0 0 0 0
8 6 1
. & 1 B 3 0
4 105 108 105 105 0 0
5 384 400 140 20 1 0
945 945 945 945 945
6 3840 4384 1800 340 30 1
10395 10395 10395 10395 10395 10395

15.7.5 Asymptotic Normality

With the recurrence relation (15.3), we can derive the asymptotic normality for X,.
Note that, X, can be expressed in terms of the sum of n independent but not
identically distributed Bernoulli random variables, i.e.,

Xn=h+1L+- -+ 1,
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where I, is a Bernoulli random variable with the distribution given by

1
P(I :1):MZI_P(ImZO)’m:LZN",n-

Therefore, we have the following asymptotic normality.

_ d
Theorem 15.1. Asn — oo, X2=E&En) % N0, 1).

N Var(Xy)
. . . _ Im—E(nm)
Proof. We consider the normalized random variable Z, ,, = oA Then
E(Zym) = 0 and anzl EZ,%,m = 1. In addition, since VarX,, = O(Inn), for

any fixed ¢ > 0, taking sufficiently large 1, we have (Inn)~'/2

that, for any fixed ¢ > 0,

< &, which implies

n
Z E(|Zn,m|2; |Zn,m| >¢g) — 0,

m=1

as n — oo. By the Lindeberg—Feller Central Limit theorem, for example, p. 116
in [3], the asymptotic normality holds.

15.8 Conclusions

There is a plethora of discrete probability problems coming from a variety of
research disciplines. We have presented a sample of these problems. We have devel-
oped a probabilistic recurrence relation methodology to solve these problems. There
are some problems remain unsolved.
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Chapter 16

On Some Inequalities

of Chernoff-Borovkov-Utev Type
for Circular Distributions

B.L.S. Prakasa Rao

Abstract We discuss some classical inequalities such as Wirtinger inequality and
weighted Wirtinger type inequality for 2 -periodic functions and study their appli-
cations for obtaining Chernoff-Borovkov—Utev type inequalities for probability
distributions with support [0, 277]. In addition we derive Chernoff type inequalities
for the wrapped normal distribution and von-Mises distribution.

16.1 Introduction

Let X be a standard normal random variable and g(-) be an absolutely continuous
function. The inequality

Var[g(X)] < E([g'(X)]?) (16.1)

was first proved by [9] and later rediscovered by [4] as a special case of a general
result for log-concave densities and was obtained independently by [5]. Analogous
inequalities for several discrete and continuous distributions are known. A matrix
variance inequality for the normal distribution was recently obtained by [10]. Matrix
variance inequalities for some multivariate distributions are derived in [13]. For a
recent overview on some stochastic inequalities and stochastic identities, see [14].
Borovkov and Utev [3] obtained a general inequality of the type (16.1). We will
discuss similar inequalities for random variables defined on the interval [0, 2r]. It
is known that the concept of mean and variance for circular random variables do
not have the same interpretation as they do for random variables taking values in
the real line. Statistical methods for analysis of directional or angular data are of
great interest. Following [6], we derive some inequalities for circular variance for
circular random variables and obtain some inequalities for standard wrapped normal
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distribution and standard von-Mises distribution. Derivation of bounds on moments,
in particular on variances, for functions of circular random variables is of interest in
directional data analysis as the exact distributions of these random variables might
be difficult to compute as in the usual case of functions of random variables tak-
ing values in the real line. The variance bounds for functions of circular random
variables derived in Sect. 16.3 deal with the general case. However it is possible
to derive better bounds for the wrapped normal distribution and von-Mises distri-
bution. These are discussed in Sects. 16.4 and 16.5 respectively. We now discuss
some classical inequalities from analysis such as Hardy’s inequalities with weights,
Wirtinger inequality and weighted Wirtinger type inequality which are also related
to the problems we are investigating.

16.1.1 Hardy’s Inequalities with Weights

Hardy’s inequality (cf. [16, p. 20]) states that if p and b satisfy the conditions 1 <
p < ooandbp < —1, then

00 p 1/p 00 » 1/p
[ / dx] < [ / 41 o) dx] (162)
0 0

T bp+1
and the constant bp_% is the best possible. Muckenhoupt [8] generalized this

inequality and proved the following result.

b * d
x/of(t)t

Theorem 16.1. Suppose . and v are Borel measures and 1 < p < oo. Then there
is a finite constant C for which

L

if and only if

D 1/p 00 1/p
du(x)} sc[/o If(x)l”dv(X)] (16.3)

1/q

_ \p r ﬂ)—t]/P
B = sup (. 0) [/0 ( @) ax (16.4)

where Z—; denotes the Radon—Nikodym derivative of the absolutely continuous com-
ponent of v with respect to the Lebesgue measure and q is such that % +1 =1

q
Furthermore, if C is the least constant for which (16.3) is true, then

B <C < p'/P1Vap (16.5)

for p>1land B =C forp = 1.
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An extensive discussion on inequalities of this type and related inequalities
known as the logarithmic Sobolev inequalities is given in [2].

The following discussion deals with inequalities for 2m-periodic absolutely
continuous functions.

16.1.2 Wirtinger Inequality

Suppose g is a 2 -periodic and absolutely continuous function with g’ € L?[0, 27)
where g’ denotes the derivative of ¢ whenever it exists. Further suppose that

2n
[ g(0)do = 0.
0

Then it is known that

2n 27
[ [g@)Pdo < [ (¢ (0)2d0
0 0

equality occurring if and only if g(6) = Acos(6 — y) for some constant A and
0 < 6y < 2x. This inequality is known as Wirtinger inequality. This can be derived
easily in the following manner.

Suppose {a,} denotes the sequence of complex Fourier coefficients of the func-
tion g. Then those of g’ are {ina, }. Applying Parseval’s identity to g’ and observing
that

1 2n
ap = —/ g(0)do = 0,
21 0
we get that

oo

1 2 oo 1 2
o [0 KO = 3 wlaal = Y lanl? =5 [0 HORA

n=—0oo n=—0oo

This result has been recently extended by [15].

16.1.3 Weighted Wirtinger Type Inequality

Let C(a, b) > 0 denote the best constant in the weighted Wirtinger type inequality

2w 2w
/0 [5(0)Pa(6)dd < C(a.b) /0 £/ (0)12b(6)d6 (16.6)

where g is a 2z -periodic function and

[mgwmwmezu
0
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Let B(L) be the class of functions a(-) € L°°([0,2m]) such that a(-) is 2x-
periodic and
inf a(@)=1and sup a(f)=L.
0<6=<2m 0<0<27
Suppose a(0) = [y(0)]? and b(0) = [y(0)]? for some y(0) € B(M),M > 1
and p,q € R such that p + g > 0. Then the weighted Wirtinger type inequality
(16.2) holds with

2 _ )
C(a,b) < |:% 0 [y ()] q)/2d9:|

2 arctan(M —(P+a)/4)
from [15]. Suppose p = 0 = ¢ and a(f) = b(A) = 1. Then it follows that

C(1,1) < 1 leading to the classical Wirtinger inequality. Suppose that b(0) = a(0)
and p = g = 1. Then it follows that

2
C(a,a) < JIT—6[arctan(L_1/2)]2

proved in [11].

16.2 Borovkov-Utev Inequality

Suppose V¥ is a random variable with a distribution function
V(@) =aVi(6)+ (1 —a)l2(0),0 <0 <2n

where 0 < o < 1 and further suppose that V; has the density function v{(6). In
addition, suppose that, for some 6y in [0, 27] and ¢ > 0,

21
[ (0 — 60)V(d0) < evi (@), by < ¢ < 27 167
[
and ’
[ (B0 — O)V(d8) < evi(@), 0 < b < b. (16.8)
0

Theorem 16.2. Suppose the conditions (16.7) and (16.8) hold. Then, for any abso-
lutely continuous function g on [0, 2],

Elg(¥) - g0 = ~E(g'W)P). (16.9)

The inequality (16.9) follows as a special case of Theorem I in [3]. We give the
proof here for completeness.
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Proof. Observe that

2 0 2
E([g(y) — g(00)]*) = [0 [ [0 g'(¢)d¢} V(d9) (16.10)

2

-/ 2 [ / 0 g’(¢)d¢} V(d6)

2

+ [O ” [ [0 ” g'(¢)d¢} V(do)

2 2]
< / [(9—%) / (g’(qs))zdﬂ V()
6o 6o
8o 8o
4 /0 |:(90 —0) /9 (g/(qs))zdﬂ V(do)
2w 2w
_ [9 [(g/(m)z [ <9—90>V(d9>] d¢
6o ¢
+ /0 [(g/(m)z /0 (o —9)V<d9)} dp
21 90
<o [ @@Pn@as+c [0 (¢ (#)°71(9)dg

2w
=<, (&' (@)*vi(p)de

=

E([g'()P).

RIS

The above inequality also holds for distribution functions on R with support not
necessarily compact under conditions similar to those in (16.7) and (16.8) (cf. [3]).
As a particular case of Theorem 16.1, the following result can be obtained. O

Theorem 16.3. Suppose ¥ is a random variable with density function v(-) on
[0,27] and there exists 8y in [0, 2] and ¢ > O such that

2w

(6 — Bo)v(0)dB < cv(¢), B < ¢ <27 (16.11)

and ;
/ (B0 — 0)v(0)dO < cv(¢h),0 < ¢ < . (16.12)
0

Then, for any absolutely continuous function g(-) on [0, 27],

Elg(¥) — g(60)]* < c E([g'(¥)]). (16.13)
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The following result follows from inequalities derived in [3] following the
literature on differential equations.

Theorem 16.4. Suppose  is a random variable with the density function v(-) on
[0,27] and there exists ¢ > O such that either

2r
g(0) = 0,/ v(p)dp <cv(0),0 <6 <2xn (16.14)
[%
or ;
gn) = 0,/ v(g)de < cv(0),0 <0 <2xm (16.15)
0

where g(+) is an absolutely continuous function on [0, 2x]. Then

E([g()]?) < 4c*E([g (W)P). (16.16)

The following two theorems give bounds on the second moments of absolutely
continuous functions of random variables defined on the real line.

Theorem 16.5. Suppose X is a random variable with the distribution function F
and the density function f > 0 on R and g is an absolutely continuous function
with E([g'(X)]?) < oo. Further suppose that

Jim g?()F(x) = lim _g*(x)F(x) = 0.

Then there exists a positive constant Cg depending on the function g and the
distribution function F such that

(E[g2(X)])? < CoE([g' (X))

Proof. Applying integration by parts, we get that

/ g2 (x) f(x)dx = [¢%(x) F(x)]% —2/ g)g () F(x)dx  (16.17)

= _2/_ g(x)g' (x)F(x)dx
-2 SO 505 o) VT dx.
e S

Applying the Cauchy—Schwartz inequality to the integral on the right side, we get
that

00 2 0o ,2 2 00
2 [T E0F ,
[ ewsemar] < [T DT roas [ wirema

equality occurring if and only if
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g(x) F(x)

o and g'(x)
are linearly related. In other words
(E[g2(X))? < CLE([g' (X)) (16.18)
where X F(X)
e =45 (S5
O

Remark. The inequality (16.18) is not of major interest as the constant C depends
on the function g(-).

Theorem 16.6. Suppose X is a random variable with the density function f > 0
on R and g is a continuously differentiable function with E([g(X)]2) < oco. Further
suppose that f is continuously differentiable such that

| llim f(x)gx) =0.

Then
(E[g'(X)])? < E[g>(X)II(f)

where 1(f) is the Fisher information.

Proof. Letu = \/7 and i = g. Note that 4 and / are continuously differentiable
functions such that
lim w?(x)h(x) = 0.

|x]—>o00

Benson [1] proved that (cf. [7])
oo
/ ([ () + (' (x) + h?(x))u?(x))dx = 0.
—00
Replacing i by Ak and applying the above inequality, we get that
oo o0 oo
/12/ h?(x)u?(x)dx + k/ I (x)u?(x)dx + / [ (x)]?dx >0
—00 —0o0 —00

for all A € R. Hence

) 2 00 00
U h'(x)uz(x)dx} 54[/ hz(x)uz(x)dx] U (u’(x))zdx].

Then it follows that
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00 2 00 00 ’ 2
[ ermax] <[ [~ s [ [ (Z3) f(x)dx}

or equivalently

E[g'(X))? < E[g*(X)(f)

where I( f) is the Fisher information. O

Remark. This inequality is the Cramer—Rao inequality. Other inequalities of these
types are derived in [12].

16.3 Circular Random Variables

Let Z be a random variable with values on the unit circle. We identify Z with a
random variable ¥ such that Z = eV, 0 < ¥ < 2m. For convenience in notation,
we denote the typical values taken by ¥ by 8. Let V' be the distribution function
of ¥. Then V(0) = 0 and V(27r) = 1. We extend the function V to R by the
equation V(6 + 2m) — V(6) = 1. Suppose V is absolutely continuous with the
probability density function v(-). Then:

(a) v(0) >0,—c0 < 6 < 00
(b) v(B 4+ 27) =v(0),—00 < 8 < 00
© [T v(@)do =1
(cf. [6]). Suppose there exists y such that
2w
(0 — 6p)v(0)do = 0. (16.19)
0

Then the conditions (16.11) and (16.12) hold provided
2w
/ (0 — 0p)v(0)dO < cv(¢),0 < ¢ <2m. (16.20)
¢

This can be seen by checking that

2w

¢
/ (6o — 0)v(0)do = (6 — 6p)v(0)dO (16.21)
0 ¢

for 0 < ¢ < 2m. Hence for any absolutely continuous function g(-) on [0, 27],

Elg(¥) — g(00))* < cE(Ig' ()] (16.22)

whenever (16.20) holds.
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Suppose ¥ is a circular random variable as discussed earlier with density func-
tion v(-). Then u is called the mean direction of ¥ if

Efsin(y —p)] =0

o is called the circular variance if
02 =E[l —cos(yy —p)] = E |:25in2 (W_;H)} .
We now obtain an upper bound for

ele((*5")) 0]

where g(-) is an absolutely continuous function. We follow the technique used in
proving Theorem 16.1.

Theorem 16.7. Suppose V¥ is a circular random variable with density function v on

[0,27]. Let
60— 6,
A(0) = 2sin (%) (16.23)
for 0 < 6,60y < 2m. Further suppose that there exists ¢ > 0
2w
A(O)W(0)dO < cv(t),t > Oy (16.24)
t
and ,
/ A(O)W(0)dO < cv(t),t > 6. (16.25)
0

Let g be an absolutely continuous function. Then
_ 2 _ 2
s (o(52)) o] s (254

Proof. Note that

2
E |:g (sin (‘/’ ; 9")) - g(O):| (16.27)
1 .
5 O

(5%

(16.26)
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[ 2 (5]
[ (5] o
L [ (o2
L] oo

The proof follows from the inequalities derived in (16.27) and an application of
Fubini’s theorem. |

dzv(&)d0:|

The following inequality follows as a special case from the weighted Wirtinger
inequality stated in Sect. 16.3.

Theorem 16.8. Suppose V is a circular random variable with a bounded, 27 -
probability density function f on [0,27x]. Further suppose that inf f =L > 0
and sup [ = M < oo. Then, for any absolutely continuous function g(-) which is
2 -periodic,

E(g(W)?) = C(f£. NHEIE' W) (16.28)
whenever E[g(¥)] = 0 where
2
C(f f) < %(arctan(Ll/zM_l/z))_z. (16.29)
Proof. Let a = and b = 4. Then a = b is bounded, 27 -periodic and inf, = 1

and sup, = L . By the welghted Wirtinger inequality, it follows that

21 2
/ [5(O)Pa(®)do < (a.a) / a (8¢ (6)12d6
0 0

if g is 2m-periodic and
2r
/ g(0)a(0)dd =0
0

where
2

C(a,a) < %(arctan(Ll/zM_l/z))_z.

Rewriting these inequalities in terms of the function f, we obtain the inequality
stated in (16.28). |

Remark. Ricciardi [15] proved that equality holds in (16.28) if and only if
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A

T=f(9+¢)

for some ¢ in R where

f@) = 1 if0<6<(x/2), andw <6 < (37/2)
=M/Lif (7/2) <60 <m, and 3/2) <0 <2m.  (16.30)

Furthermore equality holds in (3.10) with @ = f(0 + ¢) if and only if g(9) =
g(0 + ¢) where

2(0) = sin[ﬂ(@ — %)]ifo <6< g (16.31)
= (M/L)""? cos [ﬂ (9 — 377{)] if% <f<m

=—sin|:\/ﬁ(9—5%)]ifn <0< 377[
= —(M/L)""?cos [ﬂ (9 — 7%)] if 3771 <6 <2m
where . = (72 /16)(arctan(M/L)™?).

It is clear that the inequality (16.28) in Theorem 16.8 can be restated in terms of
the variance of the random variable g () in the form

Var[g(¥)] < C(f. /E(g W) (16.32)

under the same conditions as stated in Theorem 16.8 without the additional condi-
tion E[g(y)] = 0.

16.4 Chernoff Type Inequality for Wrapped Normal
Distribution

A circular random variable ¥ is said to have the standard wrapped normal distribu-
tion if its probability density function is of the form

v(0) = @2m)"* > exp (—%(9 + an)z) , 0<6<2m (16.33)

k=—00

This distribution is unimodal and is symmetric about & = 0. It is possible to obtain
a Chernoff type inequality for the wrapped normal distribution from the inequality
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(16.1) for the normal distribution by restricting the inequality to 2z -periodic func-
tions. However we will obtain the inequality directly.

Let g(-) be any absolutely continuous function on [0, 27]. Then g(-) is almost
everywhere differentiable on [0, 277]. Let I (A) denote the indicator function of set A.
Note that

2

2]
(/0 g’<¢>d¢) ( 116 > ¢lg’ (¢)d¢) (16.34)

( 116> glas ) (| T dlle (P

— 6 (/02”1[9 > 1l $)] d¢)

IA

Hence
0 2 2w
E ( / g’<¢>d¢) <E [9 ( [ 6> ¢][g/<¢)]2d¢)} (16.35)
0 0
2
- /0 EOI[9 > ¢)[¢ ($)Pd
Therefore
27
El(g(6) - g(0)?] < /0 E@I0 > ¢Dlg' @)Pdp.  (1636)
Observe that
o 1 )
E@OI[0 > ¢]) = J__ > / 6 exp (—5(9 + 2k7) )d9 (16.37)
_ LTy 1 2
=75 ), k;ooOexp( 2(9+2k7[) )d@
For k > 0,

2k 427

1 1
6 exp __(9+2k7{)2) do = / (o — 2km)exp (——oz2 do (16.38)
2 p+2n 2

© 1
< [ o exp (——az) da
o+2km 2

2w
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o0
oe(52)]
=|—exp(—za
2 [¢+2k7]

1
< exp (_§(¢ + 2k7r)2) .
Hence

1 2 1
— Y 0 exp (—5(9 + 2k7r)2) do  (16.39)

EOI > ¢)) = )
k=—o00

IA

J_ Z exp (——(¢ + 2km) )

1(¢p) + — \/_ Zoexp (—5(9 + 2k7r)2) (say).
Combining the inequalities (16.36) and (16.39), we get that
2w
E[(g(6) —£(0))%] < [0 1(9)[g'(9)de (16.40)
v TP exp (—1(¢ + 2n)2) a9
Var Jo k=0 2

2m
5[0 @) (@)Pdp + E([g' (D).

16.5 Chernoff Type Inequality for von-Mises Distribution

A circular random variable ¥ is said to have the von-Mises distribution if it has a
probability density function of the form

fO, k)= explk cos(60 — u)],0 < 6 <2x (16.41)

210 (k)

where k > 0 and 0 < u < 2. Here the function /o (k) is the modified Bessel
function of the first kind and order zero, that is,

oo

lo) =Y s (/)7

r=0

The parameter p is called the mean direction and the parameter k is called the
concentration parameter. Suppose that ;1 = 0. We denote the normalizing constant
in (16.41) by C and the function f(6,0, k) by f%(6) for convenience.
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Let g be any absolutely continuous function. Then the derivative g’ exists almost
everywhere. Then

0
g(sinf) — g(0) = [ g'(sinu) cosudu (16.42)
0

for0 <6 < 2. Let

h(u) = 1 if cosu > 0, (16.43)

=—1if cosu <0
for0 < u < 2m and

p@,u) = h(u)cosuif 0 <u <86 (16.44)
=0 otherwise.

Then

0 2
[g(sin ) — g(0)]* = /0 g’ (sinu) cosu dui| (16.45)

. ¥ ¢'(sinu) 2
= [0 n) h(u) cosu du

r P2 /(o
— / g g dui|
LJO

2

h(u)

r r2m 2 s 2
[0 P(9,u)du] |:/; (g}(ls(l;)u)) P(9,u)du:|

IA

2w 2r
= / p(6,u) du] |: (g’ (sinu))? p(6, u) du]
0 0

(since h(u) is either +1 or —1)

27
= )’(9)[ | (g'(sinu))? p(6,u) du] (say).

Hence

2n

E([g(siny) —g(0)]*) < E (V(I/f)[ | (¢'(sinu))*p(y, u)du]) (16.46)

2
- [0 Ely () p(y. 0)(g' (sinu))? du,

Observe that
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27
Ely(W)p(, )] = /0 y(0)p(0.u) fr(0) db (16.47)

2w
< / Y (8)h(u) cos u fi (8) 6

u

2w
= h(u) cosu/ v(0) f1(0) db.

u

Combining the inequalities (16.45) and (16.46), we get that

27 27
E((g(siny) - gO)) < [0 [h(u)cosu{ / y(@)fkw)de} (g’(sinu))z}du.

(16.48)
Note that
2w 2m t
[ y(t) fr (t)dt = fr (@) |:[ h(u) cosu du] dt (16.49)
0 0 0
2w 2w
= / |: fk(t)dt] h(u)cosudu
0 0
2w
= [ [1 — Fr(u))h(u) cosudu
0
(where Fi(0) = /00 fr()dt,0 <0 <2m)
0
2w
d
< /0 | cosuldu
<2r -6
< 2m.
The above inequalities show that
2w
E([g(siny) — g(0)]?) < 27 / | cos u| (g’ (sin u))*du (16.50)
0
< 27 E[(g'(siny))?| cos | (fx (¥)) 1.
Note that f;(6) > Cke_k forall 0 < 6 < 2x. Hence
: 2 27Tek /(s 2
E([g(siny) — g(0)]) = C—kE[(g (sin y))”| cos ¥/|]. (16.51)

Remark. The Chernoff type inequality for the von-Mises distribution can also be
derived from the classical Wirtinger inequality by the following method. Suppose
g(+) is a 2m-periodic absolutely continuous function with g’ € L?([0,2x]) and
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further suppose that

2w
/ g(0)do = 0.
0

Note that

A

Var[g(¥)] < E[g(¥)] (16.52)
27
< /O SO fi(6)d6

A

IA

2
sup fi () [0 g(6)]2d6

IA

2w
Supfk(9)/0 [¢'(0)]%do
sup /() [?
inf fx(0) Jo

sup fx(6) L2
WE(L%’ ¥)1°)

= exp(2k) E([g'(¥)I*).

[g" (O] fic(8)dO

IA

IA

In particular,

A

Var[g(sin(y))] < exp(2k) E[(g' sin(¥)))? cos*(y)] (16.53)
exp(2k) E[(g'(sin(y)))?| cos(y)]].

IA
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Chapter 17

Revisiting Local Asymptotic Normality (LAN)
and Passing on to Local Asymptotic Mixed
Normality (LAMN) and Local Asymptotic
Quadratic (LAQ) Experiments

George G. Roussas and Debasis Bhattacharya

Abstract Let Xy,..., X, be a random sample of size n from an underlying para-
metric statistical model. Then the basic statistical problem may be stated as follows:
On the basis of a random sample, whose probability law depends on a parameter 6,
discriminate between two values 6 and 6* (6 # 6*). When the parameters are suf-
ficiently far apart, any decent statistical procedure will do the job. A problem arises
when the parameter points are close together, and yet the corresponding probabil-
ity measures are substantially or even vastly different. The present paper revolves
around ways of resolving such a problem. The concepts and methodology used are
those of contiguity, Local Asymptotic Normality (LAN), Local Asymptotic Mixed
Normality (LAMN), and Local Asymptotic Quadratic (LAQ) experiments.

17.1 Introduction, Notation, and Assumptions

A brief description of the organization of this paper is as follows. In Sect. 17.1, much
of the needed notation is introduced, and assumptions under which a set of basic
results, Theorems 17.1-17.7 and Theorem 17.8, are established. Some statistical
applications, pertaining to testing hypotheses, are given in Sect. 17.3. In Sect. 17.5,
some applications of Theorem 17.8, relative to asymptotic efficiency of estimates,
are presented, whereas the next section is devoted to various comments. General-
izations of Theorems 17.1-17.7 and Theorem 17.8 are presented in Sect. 17.7. In
the next section, the concepts of LAMN and LAQ are introduced, and then a set of
assumptions, under which LAMN obtains, is given. In Sect. 17.9 some basic results
under the LAMN framework, Theorems 17.15-17.19, are given. In Sect. 17.10, two
examples, pertaining to the LAMN case, and one example pertaining to the LAQ
case, are discussed, and the paper is concluded with Sect. 17.11, where the proofs
of Theorems 17.1-17.6 are presented.

G.G. Roussas ()
Department of Statistics, University of California, Davis, CA 95616, USA
e-mail: roussas @ucdavis.edu

M.T. Wells and A. SenGupta (eds.), Advances in Directional and Linear Statistics, 253
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Throughout the paper, all limits are taken as n — 00, unless otherwise explicitly
stated. For the sake of simplicity, it will be assumed that the underlying r.v.’s are
i.i.d.; in a subsequent section (see Sect. 17.7), generalizations already available will
be mentioned. To this effect, let Xo, X1, ..., X, be i.i.d. r.v.’s defined on the prob-
ability space (X, A, Pg), 8 € ® open C RF k> 1,let A, = o(Xo, X1,...,Xn)
be the o-field induced by the r.v.’s Xo, X1...., X, and let P, 9 = Py | A, be the
restriction of Py to A,. It is assumed that Py g and Py g+ are mutually absolutely
continuous, Py g ~ Py g+, 0,0 € © (6 # 6*), and let

q(Xo ) Pog
be a specified version of the Radon—Nikodym derivative involved. Set

0;(0,0%) = ¢(X;:0,0%) = [q(X;:6,0%)]"2, (17.1)

so that ¢; (6, 0*) is square Py g-integrable, and the likelihood and log-likelihood
functions are given, respectively, by

L(@@)_dP”

—H%w0>

An(6,0%) = log L(8, 9 ).

In the following, we restrict ourselves to 6*, which are close to 6; i.e.,

hn .
O, :9+7%W1thhn — h e Rk, (17.2)
Thus, the relevant likelihood and log-likelihood functions are
dpP
La(0,6,) = =20 A,(0,6,) = log L,(6, 6,). (17.3)
dP, g

At this point, recall the definition of quadratic mean differentiability of a random
function.

Definition 17.1. Let g(-;0), 6 € ©®, be a random function defined on the prob-
ability space (X, A, P), and let fX g%(;0) dP < oco. Then g(-; 0) is said to be
differentiable in g.m. at 6 when P is employed if

7l (g0 +h) —g(0) = 1'g(:0)] > 0inqm.[P] as (0 #)h — 0,

where g(-; 0) is the derivative in q.m. of g(-; ) at 6, a k-dimensional random vector.
Alternatively,
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! [g(:0 +Ah) —g(0)] — K g(-;0)ingm.[P]as (0 <)A — 0

>|

uniformly on bounded sets of values of 4.

Assumptions

(A1) The probability measures { Py 9; 6 € @} are mutually absolutely continuous.
(A2) (i) For each 8 € O, the random function ¢g (6, 8*) (as defined in (17.1) for
j = 0) is differentiable in q.m. with respect to 8* at & when Py is
employed.
Let ¢o(f) be the derivative in q.m. of @o(8, 6*) with respect to 8* at
(0, 0) — a k-dimensional random vector. Then

(i1) ¢@o(0) is Ap x C-measurable, where C is the o-field of Borel subsets of ©.
Let I"(0) be the covariance function defined by
r(0) = 4&lgo(0)@o(9)]. (17.4)

Then
(A3) I'(0) is positive definite for every 6 € ©.
(A4) Foreach 0 € ©:

() g(Xo;0,0%) — 1in Py g-probability, as 6* — 6.
(1) ¢(Xo;0,0%)is Ay x C-measurable.

In terms of the q.m. derivatives ¢; (), define the k x 1 random vector A,(0) by
2 n
An(8) = ﬁ;ww), (17.5)

observe that, with I"(8) defined in (17.4),
W' (0)h = 4E9[h Go(0)1>. h € R,

and set |
A(h,0) = Eh’F(Q)h. (17.6)

17.2 Some Basic Results

We are now ready to state some basic results in the first part (Sects. 17.2
through 17.7) of the paper, which are the following.
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Theorem 17.1. Let 6,, L,(0,6,), A, (0), and A(h, 0) be defined by (17.2), (17.3),
(17.5), and (17.6), respectively. Then, under assumptions (Al)—(A4),

Py,
An(6,60,) — W A, (0) =25 —A(h, 0).
Theorem 17.2. In the same setting as that of Theorem 17.1,
L[An(0) | Pnp] = N(O.I'(8)),

where I'(0) is defined in (17.4).

Theorem 17.3. In the same setting as that of Theorem 17.1,
1
L[A(8,6n) | Phol = N (—Eh'F(G)h, h'F(Q)h).

An outline of the proof of these theorems will be given in Sect. 17.11. At this
point, recall one mode of the definition of contiguity.

Definition 17.2. Forn > 0, let P, and Q, be probability measures defined on the
o-field A, in the measurable space (X, .A,). Then the sequences {P,} and {Q,}
are said to be contiguous if whenever P,(A,) — 0, A, € A, then Q,(A4,) — 0,
and vice versa.

Remark 17.1. Although contiguity can be viewed as some sort of asymptotic mutual
continuity of the pairs P, and Q,, it should be emphasized, however, that it is
possible for { P, } and {0} to be contiguous without P, ~ Q, for any n; likewise,
it is possible that P, ~ Q, for all n, but { P, } and {Q,,} are not contiguous. These
situations are illustrated by concrete examples. See, e.g., Examples 2.1 and 2.2,
Chap. 1, in [33] (also reprinted in a paperback form in 2008).

Proposition 17.1. Let {1} be a bounded sequence inR¥, and set 6 = 6+h* / \/n.
Then, under assumptions (Al)—(A4), the sequences { Py g} and { P, gx } are contigu-
ous; in particular, so are the sequences { P, g} and { P, g, }. An outline of the proof
will be given in Sect. 17.11.

On the basis of Theorems 17.1-17.3 and Proposition 17.1, we may establish the
following results.

Theorem 17.4. In the same setting as that of Theorem 17.1,

Pn,On

An(0,6,) — W A (0) — —A(h, 0).
Theorem 17.5. In the same setting as that of Theorem 17.1,

L[An(0.6n) | Pug,] = N (3h'T(0)h, K'T(O)h).
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Theorem 17.6. In the same setting as that of Theorem 17.1,
L[Aw(©0) | Pns,] = NI (O)h, T'(9)).

Remark 17.2. (i) Theorem 17.1 may be interpreted as follows: In the neighborhood
of 6, for large n, and in the sense of probability,

An(0,6,) ~ W A,(0) — A(h, ),
or by exponentiation,
L (0, 6,) ~ EFAn@®)=A0.0) (17.7)

Expression (17.7) suggests that, in the neighborhood of 8 and for large n, the
likelihood function behaves as if it were (approximately) an exponential family.
The statistical implications of this are immense. Roughly speaking, one could
use the exponential family on the right-hand side of (17.7) to set up optimal tests
about 6 or estimates of 6, and then establish the same properties in the context
of the original family of probability measures at the asymptotic level.

(i) The precise formulation of the approximation in (17.7) is the content of
Theorem 17.7 below.

(iii)) Theorems 17.2 and 17.6 do demonstrate the point made earlier that, in the
limit, neighboring parameter points may produce definitely distinct probability
measures. The same point is made by Theorems 17.3 and 17.5.

Theorem 17.7 (Rigorous formulation of the approximation indicated in rela-
tion (17.7)). In the same setting as that of Theorem 17.1, there exists a (suitably)
truncated version AY(0) of A, () such that:

EgEN 42 ® el pBa(h) 00,

Pug[A50) # Au(8)] — 0,
Pug, [A5(0) # Au(6)] — O,

and if
R p(A) = BB ® / EV4© 4P, 9. A€ A,
A
dR 7 *
so that Sk _ E" A"(e)_B"(h), h € RF ,

dPp g

then
I Pn.o, — Rup,ll — 0, (17.8)

or

n

sup {

P PV Rup|: h € B bounded C Rk} — 0. (17.9)
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17.3 Some Statistical Applications of Theorems 17.1-17.7

For the sake of illustration, consider first the case that @ is a subset of R, and con-
sider the problem of testing a couple of hypotheses; and then consider the case that
© is a subset of R¥ (k > 2) and test a simple hypothesis.

HOCR

Recall that in an exponential family, f(x;6) = C(O)ELOT®(x), x € R,
with Q () being strictly monotone, there are level-« UMP tests for each one of the
following hypotheses against the respective alternatives; namely,

H:0=0yp, A:0>0; H:0=0p A:0<086p;

H:0<60y A:60>6; H:0>6 A:60<6;
H:0<6i0r0>6,, A:0<06<60b, (91<92).

(a) For hypotheses (alternatives) for which there are level-« UMP tests in the
exponential family, we can construct level-o tests ¢, — based on A(6;),6;,
j = 0,1, 2, boundary points — which are AUMP level-« tests

(i.e., limsup[sup(Egwy, — Egpn; O € A)] <0)
among all tests w, such that
lim sup [sup (Egwy; 0 € H)] < a.

For example, for testing H : 8 < 6, against A : § > 6, at level «, the test
defined by:

1, An(go) > Cp
$n = ¢n (An(9o)) = \Vn, An(9o) =Cn
0, An(9o) < Cn,

where ¢, and y, are constants such that &g, ¢, = a, is an AUMP test of level a.
Again, recall that in an exponential family, with Q(0) being strictly monotone,
there are level-o UMPU tests for each one of the following hypotheses against
the respective alternatives; namely,
H:@l59592,A:9<910r9>92(91<92);
H29=90,A297590.

(b) For hypotheses (alternatives) for which there are level- UMPU tests in the
exponential family, we can construct tests ¢, —based on A(6;),0;, j =0,1,2,
boundary points — of asymptotic level of significance «, which are AUMP

(i.e., limsup [sup (Egwy — Egpn; O € A)] <0)
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among all tests w, of asymptotic level of significance o, which are asymptoti-
cally unbiased
(i.e., liminf[inf (Egwp; O € A)] > @).

For example, for testing H : 6 = 6 against A : 6 # 6 at level «, the test ¢,
defined by:

On = @n (An(6o))
1, A,(80) < ayor A,(6y) > by
0, a, =< An(QO) < b,

’

(an < by) with a, — —&4/2, bu — &2 (Where &, is the pth quantile of
N (0, I'(6p))) is an AUMPU test of asymptotic level of significance .

Remark 17.3. Actually, the tests referred to under (a) and (b) above are locally
AUMP or AUMPU, respectively, but they become globally so under the additional
(usually, easily fulfilled) assumption:

Assumption (AS)

Pn~ n .
An(0)) 25 oo if /u(6y —6,) — £oo, j =0,1,2.
(i)® CRF k> 1

For testing H : 0 = 0 against A : 6 # 6y at level «, simple tests ¢, (indicators
of certain convex sets in R¥) are constructed — based on A,(6y) — which enjoy
Wald-type asymptotically optimal properties. These properties are too complicated
to be described here in any detail, but their essence may be conveyed as follows:

(P1) The weighted average power over certain surfaces (which are ellipsoids) is
largest — within a class of competing tests.

(P2) The sup of the difference of the sup and the inf of the power over certain
surfaces (which are ellipsoids) — 0.
At this point, recall that the envelope power function of a class F of level-« test
functions ¢ is denoted by B(6;«) and is defined by B(0; ) = sup{B(9; ¢);
@ € F}, where B(0; ¢) is the power of ¢ at 6 € @. Then the next property is

(P3) The sup of the difference between the envelope power and the power over
certain surfaces (which are ellipsoids) — of the test ¢, — compared to the same
sup of any other competing test have a difference whose limsup is < 0.

The asymptotic optimality properties of the test ¢, are implicit in the formulation
of (P1)-(P3).

17.4 A Convolution Representation Theorem

In this section, we consider a class of estimates, so-called regular estimates, a nor-
malized version of which is assumed to converge weakly to a probability measure.
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Then this limiting probability measure assumes of a certain convolution representa-
tion.

Definition 17.3. With 9, = 0 + Ln, the estimate T}, is said to be regular if

L [\/E(Tn - en) | Pn,Gn] = ‘C(Q),
a probability measure.
Then the following result holds.

Theorem 17.8. In the same setting as that of Theorem 17.1, and for regular
estimates,

L(O) = N, I7H(0)) * L*(0),
where L*(0) is a specific probability measure (arising in the proof of the theorem).

Thus, the limiting measure is most concentrated when it is restricted to its normal
component, and is diffused otherwise.

There are many significant applications of this theorem. Presently, we restrict
ourselves to its use in the quest of asymptotically efficient estimates; this is done in
the following section.

17.5 Some Applications of Theorem 17.8 in the Quest
for Asymptotically Efficient Estimates

Asymptotic efficiency of an estimate is looked upon in two ways: The Weiss—
Wolfowitz approach based on the asymptotic concentration of probabilities over
certain classes of sets; and the classical approach based on the variance or covariance
of the asymptotic distribution of the estimates.

The search for an asymptotically efficient estimate may be divided into two parts:
First, establish a bound for the limit (lim sup/lim inf) of a certain desirable quantity,
defined for each estimate of a target class of estimates; and second, try to identify
a member of the target class for which this bound is attained. Presently, we restrict
ourselves to the first part only.

17.5.1 The Weiss—Wolfowitz Approach

In this framework, there will be four results presented.

Theorem 17.9. For ® C R, consider estimates T, such that P, g [ﬁ (T,,—(?)fx]
— Fr(x;0), ad.f, continuously in ® for each fixed x € R and continuously on R
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foreach 0 € ©. Let L7 (0) and ut (0) be the “smallest” and the “largest” medians
of the (continuous) d.f. Fr(-;0). Then, for every 6 € ©,

t +€T(9)§Tn§9+l_2+w(9)

NG Vnoo

lim P, ¢ (9 - ) =< B(6:n.12),

where
B(0;t1,t2) = @[t,0(0)] — @[t10(0)] forallt;,t, > 0,

@ is the d.f: of N(0,0%(0)), and 6*(0) = 4Eg [¢0(6)]* . (17.10)

(Recall that ¢ (0) is the q.m. derivative at 6 of the square root of the p.d.f. of the
underlying i.i.d. r.v.’s.)

Now, allow ® C Rk, and consider estimates 7, such as those in Theorem 17.9;
ie., Py [v/n(Ty —0) <x] — Fr(x:;6), ad.f, continuously in @ for each fixed
X € Rk, and also continuously in R¥ for each # € ®. Then it is clear that, for
each h € R¥, the Py [ﬁh’(Tn —0) < x] converges to a d.f., Fr(x;0,h), say,
continuously in @ for each fixed x € R¥, and also continuously in R¥ for each fixed
6 € ©. Furthermore, let £7(0, h) and ur (6, h) be the “smallest” and the “largest”
medians of the (continuous) d.f. Fr(-; 6, k). Then the following result holds.

Theorem 17.10. For every 6 € ® C R* and each h € R¥, in the notation just
introduced and the assumptions made, it holds

lim Py g [—t1 + £7(6,h) < /ah'(Ty — 0) < t2 + ur (6, h)]
< @lno ' (0.h)] — P[—107 (8. h)],

for all t,ty > 0, where @ is the d.f of the N(0,062(8,h)), and 0%(0,h) =
W T~1(6)h.
(Recall that I"~1(#) is the inverse of the covariance I"(#) defined in (17.4).)

Next, for estimates 7,,, where a properly normalized version converges weakly
to a probability measure which has zero as its median, a result similar to that in
Theorem 17.9 holds. More specifically, we have

Theorem 17.11. Let ©® C R, and let T, be an estimate such that L]/n(T, — 0)|
P, 0] = L1, a probability measure having 0 as its median. Then

, t t
limsup P, ¢ (9 - ﬁ <T, <0+ ﬁ) < B(0;11,12),

forall ty,t, > 0, and almost all 6 € O (with respect to Lebesgue measure), where
B(0;11,13) is defined in (17.10).

If one imposes median unbiasedness to the estimates considered, then a result
such as that of the preceding theorem also holds. That is, we have
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Theorem 17.12. Let ® C R, and let T, be median unbiased estimates; i.e., for
every 0 € © and all n,

1
Pn,H(Tn > 0) > 5 and Py(T,, < 0) >

N =

Then, forall ty,t > 0 and all 0 € O, one has

, t t
limsup P, ¢ (9 - ﬁ <T, <0+ ﬁ) < B(0;11,12),

where B(0;11,t,) is defined in (17.10).

Remark 17.4. In all three cases considered above, the desirable properties of the
estimates entertained are built into the definition of the classes of estimates. How-
ever, how the convolution Theorem 17.8 is entering in establishing the inequalities
is not easy to explain without introducing many technical details. It suffices only
to say that the concentration of probabilities under the measure N (0, "1 (#)) is at
least half as large as the respective concentration under the measure £(6). This is
the decisive point on which the proofs hinge.

17.5.2 The Classical Approach

In this subsection, we state two results, each one for the case that ® C R and
® C R¥. More precisely, we have

Theorem 17.13. Let ® C R, and let T,, be an estimate such that

(i) Either Py [ﬁ(Tn —-0) < x] — &7 (x;0) forallx e Randall 8 € O,
(ii) or Py [ﬁ(Tn —-0) < x] — @ (x; 0) continuously in 0 for each x € R, where
@7 (1 0) is the d.f. of the N(0,0%(0)), for some variance 67.(6).

Then one has, respectively,

) 0%(9) > 1/02(0) and liminf [n&g (T, — 9)2] > 1/02(0), for almost all 6
(with respect to Lebesgue measure), and
(i) 0%(0) = 1/02(0) and liminf [nE(T, — 0)*] = 1/0%(0), for every 6 € ©.

For the multiparameter case, there is the following generalization of parts of
Theorem 17.13. Namely, we have

Theorem 17.14. For6 € ® C Rk, let T,, be an estimate such that
Py [Vn(T, —6) < x] = &®(x;Cr(6))

continuously in O for each x € Rk, where ®® (-;Cr(0)) is the d.f. of the
N(0, Cr(0)) with Cr(0) positive definite. Then, with I'(0) = &g [(j)o(@)(/')(’)(G)],
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Cr(0) — I'"1(9) is positive semi-definite for almost all 0 (with respect to Lebesgue
measure). Also, C(0) is continuous on ®. Furthermore, if I'(0) is also continuous,
then C(9) — I'"1(0) is positive semi-definite for all 6 € ©.

There are conditions which ensure continuity of I"(0). To this effect, make the
following assumption.
Assumption (A6)
There exists § > 0 such that, for each 8 € ® and each & € R¥, there is a
neighborhood of 6, n(0, k), with the property

248

Eg+ < M(8,h)(< o0), forall 0* € n(6, h).

h' 9o (67)]
Then we have the following result.

Proposition 17.2. Under the assumptions (Al)—(A4) and (A6), I"(0) is continuous
on 6.

To Theorem 17.14, there is the following easy but useful corollary.

Corollary 17.1. Let g be a real-valued function defined on R¥, and suppose that
(0/00,)[g0)], j = 1,...,k exist and are continuous on ©. Estimate g(0) by S, =
g(Ty), where the estimate T, of 0 is as in the theorem. Then one has:

(i) LLVN]Sn—gO)] | Pug} = N(0.03(0)), forall 6 € O, provided o3 (6) > 0,
where 05(9) =g'(0)Cr(0)g(0) and

0 0 !
#(6) = (%gw),...,@g(e)) .

(i) 0§ 8) = g'(0)I~1(0)g(8) for almost all O (with respect to Lebesgue mea-
sure).
(iii) The inequality in part (ii) holds for all 6 € O if I'(0) is continuous on ©.

17.6 Some Comments

The results stated in the preceding sections depend heavily on LAN and contiguity.
These concepts have been central in much of Le Cam’s research work, were intro-
duced by him in the literature, and were given a definite form in his seminal paper
[24]. They also constitute an essential part of his book [25], and they are found in a
more readable version in the book [26].

The important convolution representation theorem, Theorem 17.8, was estab-
lished by [17]. A version of it was also arrived at independently and at the same
time by [18]. Results employing LAN and contiguity have been obtained by the first
author in a series of papers. Relevant developments in a Markovian framework is
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the content of the book [33,38]. Contiguity was taken a step further by [15], relating
it to the statistical invariance principle.

The central assumptions among those made for the derivation of the results stated
so far is the differentiability in q.m. of the square root of the p.d.f. of the underlying
r.v.’s. Such an assumption may hold even if the pointwise derivative does not exist.
Also, it singlehandedly replaces the Cramér-type conditions involving the existence
of third-order pointwise derivatives. The interested reader may refer to the papers
[27,28].

The precise formulation of the Wald-type properties referred to in Sect. 17.3(ii)
may be found in Chap. 6 of the book [33, 38], and, of course, in the original paper
[50]. Asymptotic efficiency of estimates in terms of concentration of probability,
briefly discussed in Sect. 17.5, has been studied extensively by [51-55].

17.7 Some Generalizations of Results Stated in Sects. 17.2
and 17.4

Many of the results stated in Sects. 17.2 and 17.4 have been generalized in several
settings and different directions.

Thus, Theorems 17.1-17.4 have been carried over to the case when the underly-
ing r.v.’s are independent but not identically distributed in [29]. Theorem 17.8 was
discussed in a Markovian framework in [46], whereas statistical applications of The-
orems 17.1-17.4 and Theorem 17.8 in the same setting were presented in [21-23].
Theorems 17.1-17.4 for Markovian processes were established originally in [31],
and some statistical applications were discussed in [32,34,35]. The same theorems
were carried over to the case of general discrete time-parameter stochastic pro-
cesses in [36]. Theorems 17.1-17.4 were also discussed in the context of continuous
time-parameter Markov processes, Lévy processes, and continuous time-parameter
diffusions and Gaussian processes with known covariance by [1]. Certain aspects
of the relation between contiguity, sample size, and parameter rates were addressed
by [2, 3]. Theorems 17.1-17.4 were derived, under a sampling scheme based on
stopping times, in [4] for Markov processes, and in [5] for general discrete time-
parameter stochastic processes. The same theorems were proved in the framework
of continuous time-parameter semi-Markov processes with finite state in [39]. The
same problem was revisited by [41], where Theorem 17.7 was also established.
In general discrete time-parameter stochastic processes, various aspects of Theo-
rems 17.1-17.4 and Theorem 17.8 were discussed by [40, 44] when stopping times
are involved, and by [42,43] for fixed sample size.
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17.8 The Local Asymptotic Mixed Normal and Local
Asymptotic Quadratic Experiments

It must have become apparent by now that LAN is a valuable tool in discussing
the asymptotic distribution of the log-likelihood functions, as well as other entities
closely related to it. This is the first step in drawing statistical inference about the
underlying parameter.

It so happens, however, that LAN does not obtain in some cases which are
not too far out of the way. Two such cases are the explosive autoregressive pro-
cess of first order, and the super-critical Galton—Watson branching process with
geometric offspring distribution. What is happening instead is that, under suitable
conditions, the log-likelihood function converges weakly to a Locally Asymptoti-
cally Mixed Normal (LAMN) distribution, so-termed by [19], who first arrived at
this distribution.

For the definition of this concept, let §, be a k x k positive definite matrix such
that the matrix norm of 8! tends to 0; i.e.,

18,11 — o, (17.11)
and set
0,1, (to be shortened to 6,)) = 6 + 8;1h, h € R¥, (17.12)
so that 8, € @ for all sufficiently large n. Also, set

dP, ¢
Ay (6,,0) = log —=. 17.13
n(0n, 0) 0og AP g ( )

Then, in the language of random experiments, we have

Definition 17.4. Let§,, 6, and A, (6,, 0) be givenby (17.11), (17.12), and (17.13),
respectively. Then the sequence of experiments {(X, A,, P,g); 6 € O}, n > 1,
is said to be Locally Asymptotically Mixed Normal (LAMN), if the following two
conditions are satisfied:

(i) There exists a sequence {W,(0)}, n > 1, of A,-measurable k-dimensional ran-
dom vectors, and a sequence {T,(0)},n > 1, of A,-measurable k xk symmetric
and a.s. [P, g] positive definite random matrices, such that, for every s € Rk,

1
A (6,,0) — [h’Tnl/ 2(0)W,(6) — Eh'T,, (G)h} — 0in P, g-probability.

(17.14)
(ii) There exists an a.s. [P, g] positive definite k x k symmetric random matrix
T (6), such that,

LW (0). Tu(0)]| Png s = LAW(O). T(0)]] Pyj. (17.15)
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where W(0) ~ N(0, I) and is independent of 7'(6).
By setting,

A (0) = TH2(0)W,(6) and A(6) = TV2(0)W(6). (17.16)

properties (i) and (ii) become as follows:
() Forevery h € R¥,

An(6.0) — [ Ap(8) — ' T, (9)h] — 0 in P, g-probability,  (17.17)

(ii")
L{[AR(0). T (0)]| Pno} = LLUA®), T (0)]]Pg)}- (17.18)
If conditions (i) and (ii) (or (i’) and (ii")) are satisfied, we also say that the sequence

of families of probability measures { P, g;6 € ©}, n > 1, has the LAMN property
ateach 0 € ©.

Remark 17.5. Comparing the convergencesin Theorem 17.1 and in relation (17.17),
it is seen that the constant A(h, 0) is replaced in (17.17) by the r.v. %h’Tn(G)h.
Also, comparing the convergences in Theorem 17.2 and in relation (17.12), it is
seen that the random vector A, () is replaced in (17.18) by the random vector
[A4,(0), T,,(0)], whose asymptotic distribution, under P, g, s L{{A(0), T(0)] | Pg}
rather than N (0, I"(0)). See also Remark 17.7 for some additional comments.

Remark 17.6. There exist statistical experiments (to be discussed in Sect. 17.10,
Example 17.3), defined by (17.17) and (17.18), but for which the relations stated in
(17.16) may not hold. Such experiments are called Locally Asymptotic Quadratic
(LAQ) experiments [20, 26]. The points at which LAN or LAMN do not hold
are called ‘critical points’. For LAQ experiments, it is observed that, under the
contiguity of { P, ¢} and { P, g, }, the following relation holds:

Elexp(h’ A= LW'Th)] =1 forallh, (17.19)

where A and T are as they appear in (17.18).

The assumptions made below refer to the segment of r.v’s Xi,..., X, com-
ing from a general (discrete-time parameter) stochastic process {X,}, n > 1; this
process need not even be stationary, either in the strict or in the wide sense. In
the present context, the o-fields A, are induced in X by the segment of r.v.’s
X1, Xy

Assumptions Pertaining to the LAMN Case

A set of assumptions will be listed below which imply the LAMN property, after
some additional notation is introduced. First, it is assumed that, for j > 1, a
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regular conditional probability measure of the distribution of X;, given X; | =
(X1,...,X 1), is absolutely continuous with respect to a o-finite measure y ; with
corresponding conditional p.d.f. f;(- | X;_1; 6), and the distribution of X is abso-
lutely continuous with respect to a o-finite measure p with corresponding p.d.f.
f1(-; 8). Furthermore, it is assumed that the support of all p.d.f.’s are independent
of § € ®. These assumptions imply that, for any 8, 0* € @, P, ¢ and P, g+ are
mutually absolutely continuous, P, g ~ P, g+, n > 1.
For notational convenience, set

X 1 X-1:0) = f;(0), =2 J1(X1 | Xo;0) = f1(X1:0) = f1(0).

Assumptions
(B1) With 6, = 6 4+ ;' h and f;(6) = f;(X,|X,_1:6), define &, (-; 6, h) by

Enj (0.0 = £]200) = £]720) = £}2C1Xj20:00) = £ C 1 X000).

Then assume that there exists a k-dimensional random vector &;(6), such that

— 0,

2
[ [geeon = 5w5150) | an,

k
2 Eo
j=1

(B2) Setn;(8) =§; (9)/fj1/2(9),j > 1. Then assume that
Eolnj (@) | Aj—1]=0 as[Ps]. j =1

(B3) There exists a k xk symmetric, a.s. [ Pg] positive definite random matrix 7(6),
such that

n
5,1 € [nj(0)m;(6) | Aj—1] ¢ 8," = T(6) — 0in Py-probability.
j=1
(B4) For every ¢ > 0 and every h € R¥,
- 2
Y & {|h'3;1nj(9)| I[|W's; n;6)| > 8]} 0.
j=1
where, of course, /(-) stands for the indicator function.

(B5) Either for every h € Rk, there exists k > 0, such that
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z 2
-1
supZ&; W8, 0 (0)|" <,
n>1 i=1

or
nEy [1'8; " n; (t9)|2 = O(1), uniformly in j.

(B6) Define the 1.v.’s 1, (0, h) by:

129
@y = L0y

117%0)
Then assume that, for every h € RF ,
néy {[nnj(G,h) +1]* - 1} = O(1), uniformlyin j > 1.
The main results to be stated in the next section also require a specification of

the (fixed) matrix d,, the random matrices 7,(6), and the random vectors W, (0).
Namely, 6,, T,(0), and W,,(0) are taken to be as follows:

n

Spn =Y € [n; ()0 (6)]. (17.20)
j=1
Ta(0) = 8," 3> " Eo [0y (O)n;(0) | Aj—1] ¢ 6, (17.21)
j=1
Wa(0) = T,7'2(0) | 8, D " n;(6) | - (17.22)
j=1

17.9 Some Basic Results

Here is the first result.

Theorem 17.15. For every h € R¥, let 6, be defined by (17.12), and let 8,, T (6),
and W, (0) be defined by (17.20), (17.21), and (17.22), respectively. Then, under
assumptions (BI1)—(B6), the sequence of families of probability measures { P, g; 6 €
®}, n > 1, has the LAMN property, that is,

Ay (6y,0) — [h’Tnl/z(O) W, (0) — %h/Tn (G)h] — 0in P, g-probability for every

h e RF, (17.23)

and



17 Revisiting LAN and Passing on to LAMN and LAQ Experiments 269
LW (0), Ta(O)] | Ppot = LUW.T(0)] | Po} . (17.24)
where T(0) is as in assumption (B3), W ~ N(0, I) and is independent of T (9).

Furthermore,

Theorem 17.16. In the setting of Theorem 17.15, it holds
L[An(0n.0) | Pug] = L [h’Tl/z(G)W — LW (O)h | Pg] ,
and therefore
c [h'T,}ﬂ(e)W,,(e) — LW T, (0)h | P,,,e] Ny [h’Tl/z(G)W — LY (6)h | Pg] .

The following is a contiguity result. Namely,

Proposition 17.3. In the setting of Theorem 17.15, the sequences of probability
measures { Py g} and { Py, g,}, n > 1, are contiguous.

Theorem 17.15 and Proposition 17.3 immediately yield the following result.
Theorem 17.17. In the setting of Theorem 17.15 and Proposition 17.3, it holds
A (6, 0) — [h/T,,l/Z((?)Wn ©) - 11T, (e)h] — 0 in Py, -probability.
In the following, a certain local approximation of the underlying family of prob-
ability measures is discussed. To this end, consider A, () and construct a certain
truncated version of it, call it A% (8), like so: For 0 < k, 1 oo, define W,”

(= Wk (6)) by

W = Wal (|7,2W,

< kn) = Wal (180 < ),
and take
A T2k — T2, T (| An| < kn) = Anl (|An] < kn) .
Then it is seen that
A¥(0) — An(0) — 0in P, p-probability, (17.25)

and
Chn(0) = / exp [h/AZ(G) — %h’T,,(@)h] dP,p < o0. (17.26)
X

Next, define the probability measures

Oni(A) = Con(0) / exp [ AX(0) — LW T(0)h] dPyg, A€ A,, (17.27)
A
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so that
d Qn,h

dP, e

Then the following results may be established.

= Cpn(0) exp [ Ax(0) — ' Tw(0)h]. (17.28)

Theorem 17.18. In the notation introduced above and under assumptions (B1)—
(B6):

(@) sup[||P, g 151y — Onnll: h € B, a bounded subset of R¥] — 0.
(b) sup[|Cy (8) —1]; |h| < b] — O foreveryb > 0.
(c) ||Pn,0+b’;‘h,, — On.h, || = O for every bounded sequence {h,}, n > 1, in Rk,

It is in the sense of Theorem 17.18 (parts (a) or (c¢)) that the probability measures
P, gys-1p are approximated locally (and in the sup norm mode) by the probability
measures O j defined in (17.27). Although in the construction of the measures
On.p it was A} (6) rather than A,(6) which was used, this would be irrelevant
asymptotically, because of (17.25). The usage of A (6) was dictated in order to

ensure finiteness of the norming constant defined in (17.26).

Remark 17.7. Unlike Theorem 17.7 (see relations (17.8) and (17.9)), the approx-
imation provided here in Theorem 17.18 is not by a standard exponential family.
Instead, the approximating family is a curved exponential family, so termed by [14].
Roughly speaking, an exponential family is curved when the dimensionality of the
sufficient statistic for 6 is larger than the dimensionality of 6. For example, the nor-
mal family N (6, 62), 6 € R, is a curved exponential family. For more information
the interested reader is referred to the book [12], and in particular Sect. 16.9.

In Theorem 17.16, the limiting distribution of the log-likelihood is a curved
exponential family. However, independence of W and T'(f) and the fact that
W ~ N(0, I1) turns the limiting distribution into a normal distribution, conditioned
upon T'(6) = t. This fact is exploited for making statistical inference in the limit,
and then transposing it to the original family; optimal properties of statistical pro-
cedures will be of the asymptotic variety. Relevant references in this respect are
[6,7,13,16,47-49]. The proofs of Theorems 17.15-17.18, when stopping times are
involved, can be found in [8—11]. A thorough discussion of this sort of asymptotic
results with applications to time series models can be found in [20].

A convolution representation result similar to that in Theorem 17.8 is established
here, too. More precisely, one has

Theorem 17.19. Suppose assumptions (BI1)—(B6) hold. Also, let {V,}, n > 1, be
a sequence of k-dimensional random vectors of regular estimates of 6; that is, for
every h € R, there is a k-dimensional random vector V(0), such that

L0 (Ve = 60). Ta(O)] | Pug,} = LUAV(O). T(O)] | Po} = L(6). say. (17.29)

Let L1 gy be a regular version of the conditional distribution of V(0), given T (0).
Then



17 Revisiting LAN and Passing on to LAMN and LAQ Experiments 271

LV(0)|T(0) = LT(g) = L1 * L2 a.s. [Pe], (17.30)

where L1 = N(0, T~1(0)) and L, is the conditional distribution (under Py) of
V(0)—T~Y2(0)Z(0), given T(0), and Z(0) follows the N(0, Iy) distribution and
is independent of T ().

It is to be noted that the convolution representation here refers to the conditional
distribution of V(6), given T'(0), unlike the case in Theorem 17.8. This is the way
one may circumvent the curvature of the limiting family in the present context. A
version of Theorem 17.19, when stopping times are involved, can be found in [45].

17.10 Examples Pertaining to the LAMN and LAQ Cases

Here are three examples whose discussion falls into the LAMN and LAQ frame-
works.

Example 17.1 (Explosive autoregressive process of first order). Such a process
consists of r.v.’s X ;, j > 0, generated as follows:

Xj=9Xj_1+8j, Xo =0, |9|>1,

where the ¢ ’s are independent r.v.’s distributed as N (0, 1). The X ;’s form a Markov
process with transition p.d.f. that of N(6x,_1, 1), so that

1
N2

In [8], it has been checked that assumptions (B1)—(B6) hold, so that the underlying
family of probability measures is LAMN (see also [16, p. 110], and [49, pp. 135-
136]).

The key quantities here are:

fi(xjlxj-1:0) = f(xjlx;-1:0) = exp [—%(Xj - 9xj—1)2} :

o2 (62 — 1)h
St = TR sothat9n=9+9—n,
£y = L1O)
e
where
. 0 Xi—1(x; —0x;_ 1
fi0) = @f(xﬂxj—l;@) = %=l i/ﬂ j=1) exp |:_§(Xj —Oxj_l)z} ,

so that
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£0)  f50)

n;j(0) = fjl/z(e) =70 =xj_1(x; —0x;_1).
It follows that
ro = Co L Af W) = Zim il
(5o x2y)
An(0) = @ _I)ZX, 1€

Furthermore, it is seen that the log-likelihood is given by

(9 —Dh Yo Xj- I(Xj —0Xj-1)  (0°=1)*h? 2,
Xi_,+op, ,(1).
on (Z/=1 2 )1/2 202n ; Jj—1 .60

From results found in [6, pp. 164-165] and [16, p. 110], it is seen that

LITu(0)|Pag] = LIT(0)Po] = 17
L{AO), Ta(O)]| Pa g} = LUT > (O)W. T (6)]| P},

where W ~ N(0, 1), T'/2(9) ~ N(0,1), and T'/2(#) and W are independent (all
under Py).

Example 17.2 (Super-critical Galton—Watson branching process with geometric
offspring distribution). This example has been discussed in [6, p. 163]; [13, pp. 853—
8547; [8]; and [49, pp. 133—135]. In the present case,

1 Xj—=Xj—1 1 Xj—1
_fj(xj-|xj_1;9)=f(xj|xj_1;9)=(1—5) (5) y 0> 1.

Here, the geometric offspring distribution is given by
PXi=j)=0"'a-0"YH "1 j=1.2.. 6>1I,

so that
EgX; = 0 and Varg(X,) = 0(0 —1).

The key quantities here are (see [13, pp. 853—-854] and [49, pp. 133-135]):

5_1 _ 91/2(9 - 1)

0126 — Dh
n gn/2

, sothat 6, = 0 + on/2 )
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/i (0) 9 8 o
£;(0) = fjlj/2(9)7 where ﬁlog S(xjlxj-1:0) = log £i(6) = /9(9—_;)1’
and f;(6) = f3(6) x 55 10 f7(6), so thatn; (6) = X;— 60X
Jj J gJi nj 50— 1)

It follows that

0

1™ S (X, —0X,-
ONLES S P v 1 7Rt 721
j=1

66 —1) Gy Xj—]

and "
Zj:l (Xj - 9Xj—1)

An(e) = 9(}1—1)/2

Furthermore, the log-likelihood function is given by

h (6 —1)2h?
An(0,0) = o 1)/ZZ‘(XJ 0X ;- 1)—2—2)(, 1+ 0p, 4(1).

j=1
It can be seen (from the references just cited) that
L[Tx(0)| Pnp] = LIT ()],
L{ARO). TaO)]| Pug} = LUT 2 (O)W. T(6)]] Py},
LIAn(O)|Pap] = LITVO)W | Py,
where 7'(0) is an exponentially distributed random variable (under Pg) with unit
mean, W ~ N(0, I), and T'/2(#) and W are independent.

Example 17.3. Let us consider the process described in Example 17.1 again, where
6 = 1is the true value of the parameter 6 (a unit root autoregressive process of order
one). We use (17.20) to find §,, as indicated below, and, without loss of generality,
take 8, = n,sothat6, = 6 + %

n
> B nn—1) 2
Soxz, | ="l
j=1
We find that the log-likelihood function is given by
An(6n,0) = ZX] 1€/~ 5 ZX] L +op, ,(1).

It can be seen that the distribution of A (6,, 0) tends weakly to the limit distribution
of Ay, where
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1 h2 1 1 h2 1
A,,:[ W,th——/ Wtzdtz—(le—l)——/ W2 dt,
2 2 2
0 0 0

and W; is a Weiner process or standard Brownian motion [16,30,48]. Here

1 & 1 1
B (@) = Y Xy = /0 W, W, = SOV = 1),
i=1

where W,2 is a N(0,¢t) r.v. for fixed t,
1 ¢ 2 ! 2
T6) = 5 X7, :>/0 W2 dr,
j=1
and
1 1
L[An(0), Ta(O)] | Pnot = LI(A,T) | Pyl = (/ W, dW;, / w2 df)~
0 0

It can also be seen that

L@, = 1) | Pag) = L[3W2=1) [ [y w2 ar | Py

where ;
0= Zj:l XjXj-1
==l - -
2j=1 X7
and
Ly X (X;—Xjm1) A3 X e A (O
n(/é =1 j=147-114] =) w2j=14j-18j  Au(0)
a—1) =

Yo X7 w i Xi Ta®)

17.11 Outline of Proofs of Some of the Basic Results

From Sect. 17.1, recall that ¢ ; (6, 6*) = [q(X;:6,6%)]'/2. Keep 6 fixed, replace 6*
by 6, = 6 + hy,//n with h,, — h € R¥, and set

onj (0) = 0} (0.60) = [q(X;:0.60)]'.
Also, recall that ¢o(6) is the q.m. derivative of ¢o(6, 8*) with respect to 6* at 0

when Py is employed. In this notation and always under assumptions (A1)—(A4),
we have
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Lemma 17.1.

n

Z[%j (0) — 117 = &E[h' ¢o(0)]? in Pg-probability.
=0

Proof. See Lemma 5.1 in [33,38]. O
Lemma 17.2. With A, (6, 60,) = A, (0) defined by (17.3), we have

n n
An(0)—2 Z[gonj @) -1+ Z[(pnj (0) — 1]> = 0 in Py-probability.
Jj=0 j=0

Proof. See Lemma 5.3 in [33,38]. O

From Lemmas 17.1 and 17.2 we have, by subtraction
n
Ay (0)—2 Z[(pnj (0) — 1] = —Eg[h' ¢o(0)]* in Pg-probability. (17.31)
j=0

Now, in the alternative definition of q.m. differentiability (see Definition 17.1),
replace 6* by 6 + Jiﬁ to obtain

h q.m.
0,0+ —]—-1 Wei(0
\/E|:(pj(’ +\/’7) :|E; (pj( )»

since ¢;(6,0) = 1; or

7 [0nj (6) — 1] (%"’; 15 (6). (17.32)

4

However,
Lemma 17.3. /n[py;(6) — 1] — 2h'¢; () in the first mean [Py).
Proof. See Lemma 5.4 in [33,38]. O

Butif U, ﬂi U, then
|EUn = EVU| = |EWUn = U)| < E|Un = U| < EV2|U, —U> > 0
(see, e.g., Corollary to Theorem 2, Chapter 6, in [37]). Accordingly,
€5 [V (¢3;(0) — 1)] — E5[2h'¢; (6]

However, Sg(p,%j (6) = 1, so that E[h'¢;(6)] = 0; Thus, we have the following
lemma.
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Lemma 17.4. Foreach h € R¥, & [¢;(6)] =0.

Next, omit 6 in the expressions for ¢,;(0) and ¢; (6), and set

1 .
Y =(gnj—1)— ﬁh ©; — (Egpnj — 1),

so that £gY; = 0, and apply Tchebichev’s inequality to get the result.

IA

n
Po[ D Y| >e
j=0 i

1 - n+1 1.
8—2092 Y= 8—2092 |:((/)n0 -)- —h,%]
j=0

1
Eo |/ (gno — 1) = W go|* —> 0,

IA

so that

1 n
Z Y; = Z Onj — 1) — 7 > W¢; —(n+1)(Egpno — 1) — 0in Py-probability.

j=0 j=0

(17.33)
Combining (17.31) and (17.33), we get

Ay — «/ﬁ Z Rg;—2m+1)(Eopno—1) —> —Eo (W (po) in Pg-probability.
Jj=0

(17.34)
From the identity

Oro—1=(ono— 1> +2(pno— 1),
we obtain, by taking expectations,

0==Eg(prg—1) =Eo (9no — 1> +2(Eopno — 1), or
Eo [/ (@no — D]* + 21 (Egpno — 1) = 0. (17.35)
Since &y [ﬁ((p,,o - 1)]2 — &y (h/gbo)z, relation (17.35) yields
21 (Egpno — 1) > —Eq (Hg0)”

or .

2(n 4+ 1) (Eopno — 1) — —Eg (W' g0)” . (17.36)

Relations (17.34) and (17.36) yield then

Ap — «/_ Z Woj — —2E (W (/)0) in Py-probability. (17.37)
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Since, by relations (17.6) and (17.4),
1 1
—A(h.6) = — W T(O)h = — 2 x 48 [60(6)]” = ~285 [1'60(0)].
and
2 n
T 9O = 4O
by (17.5), then relation (17.37) establishes Theorem 17.1; i.e.,
Proof of Theorem 17.1.

An(0) —h' Ay(0) —> —A(h. 6) in P, g-probability.

We proceed now with the proof of Theorem 17.2.

Proof of Theorem 17.2. By dropping 6 for convenience, it suffices to show that, for
all h € R¥,
L (W Ap | Pyg) = N, h'Th).

From (17.5),
2 n
’ _ >
Jj=0
withther.v’s W'¢;, j =0,1,...,n,beingiid., E (h'¢o) = 0 and og (W' ¢o) < o0.
Then the CLT applies and gives

% Z Wej = N (O, &y (h’(/')o)z) under Py,
j=0

or
LK An(8) | Ppg] = N(O, K'T(9)h) forall h € R,
so that
L[An(0) | Pl = N(O, I'(0)).

Proof of Theorem 17.3. It is an immediate consequence of Theorems 17.1 and 17.2.
O
At this point, the contiguity result stated in Proposition 17.1 is established.

Proof of Proposition 17.1. By Theorem 17.3,

02 2
L(An| Ppg)= N (—7, o )
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where 02 = h'I"(9)h, and

A o’ 2 1 A o\’ 2
E*dN|(——, o =/ E* ex —()L—}——) 20 di
[l; ( 2 ) R V270 P 2 /

— 1 o\, 2 —
_/Rmaexpg—(k—T) /20§ dA =1.

Then statement (S3) on page 11 in [33, 38] implies statement (S;), which is the
required contiguity. O
Proof of Theorem 17.4. Contiguity of { P, ¢} and { P, g, } implies that, for any A, -
measurable r.v.’s T, the convergence T, — 0 in P, g-probability implies 7, — 0
in P, g, -probability, and vice versa. This is so by Proposition 2.1, page 8, in [33,38].
Applying this for A, (0) = A, (6, 8,), we obtain the desired result. |
Proof of Theorem 17.5. 1t follows by Theorem 17.3, the contiguity of the sequences
{Pn,0} and { P, g,}, and Corollary 7.2, page 35 in [33,38]. O
Proof of Theorem 17.6. It follows by Theorem 7.2, page 38 in [33, 38], by way of
Theorems 17.1, 17.2, and Proposition 17.1. O

The remaining main results are considerably more complicated, and any attempt
to provide even an outline of their proofs would take us too far afield. So, this section
is concluded here. Interested readers are referred to the different papers written by
the authors for the omitted proofs of results stated herein.
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Chapter 18
Long Range Dependence in Third Order
for Non-Gaussian Time Series

Gyorgy Terdik

Abstract The object of this paper is to define the long-range dependence (LRD) for
a Non-Gaussian time series in third order and to investigate the third order properties
of some well known long-range dependent series. We define the third order LRD in
terms of the third order cumulants and of the bispectrum. The definition of the third
order LRD is given in polar coordinates.

18.1 Introduction

Higher order spectra and in particular bispectra are very efficient tools for time series
analysis, the development of the rigorous theory has been started in the sixties [5, 8],
the applications include testing linearity and Gaussianity [14,31] parameter estima-
tions [6, 19] and so on with a huge literature. At the same time many papers have
been devoted to the research on the long memory time series. The memory of a time
series is defined usually by the order of decay of the covariance function at infin-
ity. The classical stationary ARMA models have short memory with exponentially
decreasing covariance function. The long memory means that the decay is hyper-
bolic. The asymptotic distribution of the sums of the long memory processes is well
studied due to [9, 10,23-27,32]. The dependence structure of a time series charac-
terized by the covariances only for a Gaussian series, in this case being uncorrelated
and independent is equivalent, hence long memory and long-range dependence are
synonymous concepts in time series analysis. If the time series is not Gaussian
all the higher order cumulants are necessary for the description of the dependence
structure. The independence, for instance, implies and implied by that each higher
order cumulant is zero, except all variables in the cumulant are the same. Although
Non-Gaussianity and long-range dependence have been observed in many areas, in
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network traffic [22,29,37] in asset returns and exchange rate data [11], musics [7],
etc., there are no results neither on the higher order statistics nor on higher order
spectra of non-Gaussian and long-range dependent time series.

The object of this paper is defining the long-range dependence (LRD) of a time
series in third order and investigating the third order properties of some well known
long-range dependent non-Gaussian series. We define the third order LRD in terms
of the third order cumulants and of the bispectrum. Both the third order cumulants
and the bispectrum are symmetric, therefore we consider their values on the princi-
pal domains only. The definition of the third order LRD is given in polar coordinates,
since the origin and the x-axis have some particular importance. Besides the pri-
mary singularity of the bispectrum at the origin, the singularity on the whole x-axis
is allowed. Similarly, not only the radial decay of the third order cumulants at infin-
ity is considered but its behavior when it is approaching the x-axis on the ‘circle
with infinite radius’ as well. The marginal bispectrum and the third order cumu-
lants on the x-axis couple these properties of the bispectrum and the third order
cumulants. In Sect. 18.3 we consider five basic non-Gaussian models; the fraction-
ally integrated noise, the linear fractional noise, H,-process, Rosenblatt process and
ALISDLG process serve as prototypes for the third order LRD. In the last section
we summarize the results and put some conjectures for further subjects of investi-
gations. The proofs are technical and omitted, interested readers may consult with
[36] and the author.

18.2 Long-Range Dependence

It is a well known that for an i.i.d. series X1, X5,... X, (with mean zero and
variance Var (Xy) = 0)2() the variance of the sum of Xj’s changes linearly with n,

ie.,
n
Var (Z Xk) =no%.
k=1

Asymptotically the above result holds even when Xy is a stationary time series. Let
X/ be a zero mean stationary time series given in spectral representation

12
X, = [ / elZn[a)MX (da)),
—-1/2

where E | My (da))|2 = S5 (w) dw, and S, (w) is its spectral density function. The
variance of the sum ZZ;(I) X is calculated directly from the spectral representation,
we have
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n—1 1/2 ,i2nne _
Var Z X[ = Var / WMX (da))
(=0 —1/2
1/2 2
= / Ss (w)dw.
—1/2
: : 14 i2nnw i2nw 2
Now, as far as S5 (0) is finite the Fejér kernel |(e — 1) / (e — 1)| guar-
anties the limit
1 1/2
lim — [
n—-oo n —-1/2

hence for large n we obtain

n—1
Var (Z Xe) ~nS;, (0).

=0

ezznnw _

ei2row _ |

2
S (w)dw = S5 (0),

elZnnw _

ei27rw —1

If this is true for a particular time series then it is expected that the estimated vari-
ances of the aggregated series for different n will show this approximation. Namely
estimating and plotting the logarithm of these variances against log (n) we obtain a
linear function with slope 1. Several phenomena have been reported in the literature,
see [2,28], where the slope of log (n) is different from one, usually it is greater than
1, i.e., for large n

n
log Var (Z Xk) ~ 2H log (n) + const.,
k=1

where H € (1/2,1). Half-slope H is called the Hurst exponent, the name goes
back to [17]. From now on we shall use a shifted version h = H — 1/2 of H,
since it is frequently used in time series analysis. When the slope is larger than
one, the spectrum has singularity at zero, which is equivalent to the divergence of
the covariance series Z?:—oo Cov (X¢, X¢+x)- Therefore the decay of the covari-
ances Cov (X, X¢1%) is slower, hence the dependence of the values X; and X,z
is stronger then in a usual case when the S> (0) is finite. A stationary time series X,
£ =0,+1,4£2... £ n will be called long-range dependent (LRD) if its spectrum
S> (w) behaves like |a)|_2h at zero, more precisely

S> (w)

lim ————— =qj, (18.1)
=0 o L (1/ |wl)

where & € (0,1/2) and L (-) is a slowly varying function at infinity. This definition
of long-range dependence can be stated in terms of autocorrelation function as well,
since, as far as L (-) is quasi-monoton slowly varying, (18.1) is equivalent to:
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Cov (Xe4k, Xe) _

ki)rr;o |k|2h_l Lo de, he(0,1/2). (18.2)
Note here the connection between these two constants g and g, namely the ratio
as/ae = 27272 (2h) cos (7rh) is constant. From now on this ratio will be
denoted by y,j. In other words, the autocorrelation function decays hyperbolically.
In fact although the spectrum is in L its Fourier coefficients Cov (X, X% ) are not
in L; any more. The equivalence of (18.1) and (18.2) is studied in the theory of
regular varying functions in details, we quote here the results of two Theorems,
4.3.2 and 4.10.1 of [3]. Let L (-) be quasi-monotone slowly varying function, then
forh € (0,1/2)

(o)
D L(k) [kt ek = 21 2hg=2h (21 cos () || L (1/ o))
k=—00
(18.3)
when @ — 0. The left hand side converges to a function, S» (@) say, such that it
fulfils (18.1). The Tauberian conversion of this Abelian result is also true, namely
if the Fourier coefficients of the right hand side, Cov (Xy, Xj) say, decreasing then
it fulfils (18.2). These results of calculus covers totally the problem of LRD for
Gaussian time series. It should be emphasized that either we are given the spectrum
around zero
Sx(w) =~ coyan lw| L (1) o)), w—0,

where ¢ is some (positive) constant which might depend on /4, or the covariances
at large k
Cov(Xpik. Xo) ~ co k" VL (k|), k — oo,

they defines each other including the exponents, constants and slowly varying
functions.

If the time series is not Gaussian then the higher order cumulants and spectra
provide some additional information on the structure of dependence.

18.2.1 Bispectrum and Cumulants

Let the process Xy be centered and stationary in third order, then its third order
cumulants are

Cum (X6+k1 s Xttks s Xe) =EX¢yr, Xy, Xe
= C3 (k1:2), ki,ky =0,£1,£2,...

(notation: k1.2 = (ky1,k2)). The third order cumulants are called bicovariance as
well. An easy consequence of this definition is that the following properties are
fulfilled
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Cs (k1:2) = C3 (ka, k1)
= C3(—ki,ky —k1).

These equations provide the symmetry of the third order cumulants, the plain is
divided into six equivalent parts, each of them is sufficient for the determining of
the third order cumulants on the whole plane. One of these parts, called principal
domain for the third order cumulants, is 0 < k, < kj, i.e., the lower half of the right
upper quarter of the lattice with integer coordinates in the plain.

The bispectrum S3 is a complex valued function of two variables with Fourier
coefficients C3 (k1:2), i.e.,

1/2
Cs (ki) = /] ei27r(w1k1+w2k2)S3 (1:2) dwr:s.

—1/2

In case the series C3 (k1, k») is in L; then the bispectrum has the series expansion

oo oo

Ss(@ia)= Y, Y Cs(kip)e Prekiteka) (18.4)

k] =—00 kz =—00

While the spectrum is real and nonnegative the bispectrum is generally complex
valued and since C3 (ky:») is real, we have S3 (w1:2) = S3(—wi:2), (notation:
w1:2 = (w1, w3)). The bispectrum S3 is periodic

S3(w12) = S3 (w1 +my, w2 +mz), my,my=%1,£2,...,
and it has the following symmetry

S3 (w1:2) = S3 (w2, w1)
= 83 (w1, w3).

where w3 = —w; — w;. These symmetries imply 12 equivalent domains for the
bispectrum, see Fig. 18.1, the principal domain traditionally is the triangle A with
vertices (0,0), (1/2,0), and (1/3,1/3) (A denoted by 77 in Fig. 18.1) [15,35]. The
Fig. 18.1 shows the symmetry lines going through on the origin for the bispectrum
of a continuous time process as well.

The cumulants on the x-axis will play some particular roll later on, they are the
Fourier coefficients of the marginal bispectrum M3, i.e.,

1/2
Cum (Xg. Xo. Xo) = [ M @) do
—1/2

The marginal bispectrum is calculated directly from the bispectrum
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Twelve triangles
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E 213 <12 13 0 13 12 23 1

Fig. 18.1 12 domains of bispectrum

1/2
Ms (w) = /1/2 S (@, A) dA.

The following formula is useful for the calculation of the cumulant C3 (£1:3) of
a stationary process. When the bispectrum is given we have

Cz (Ly0) = / . eizn(€1w1+€zwz)g3 (w1:2) dwi:2,
[0,1]

since the bispectrum is symmetric, see above we split the integral into 12 parts, each
corresponds to a domain A,,, m = 1 : 12, with boundary according to the symmetry
lines, see Fig. 18.1. For instance A; denotes the lower half of the upper quarter of
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the plane, it corresponds to the triangle 77 in the Fig. 18.1. Now, the transformation
from A,, into A does not change the bispectrum (conjugates it at most) it changes
the exponent only. Let us consider the integral

/ el2mtiertbam) gu (0)).5) dwya,
As

and transform As into Ay, i.e., change the variables w; = —A1 — A3, and w, = A1,
we obtain

[ e!2mlrerthm) g1 (p1,) dwry = / e 2rlmtilti=tid2) 53 (h1.5) d A2
As A
=c3(lr =41, —41),

where
s (61;2) _ / ei27r(51w1 +52w2)S3 (w1:2) dwy.
AN

After transforming all domains into the A; we obtain the following expression.

Lemma 18.1. The continuous time cumulant function can be calculated in terms of
integral c3, namely

C3(£y,£2) = 4Re (Sym ez (@1, €2) +c3 (b — L1, —41) +c3 (L1, 2 — 51)]>,

L2

where symy  denotes the sum according to all possible permutations of {1:
divided by the number of the terms.

18.2.2 Long-Range Dependence in Third Order

The relations (18.1) and (18.2) concern on the behavior of the covariances at infinity
and the spectrum at zero. Similar results in 2D are available for isotropic case only,
see [30, Ch. VII, Theorem 2.17].

Lemma 18.2. Suppose that the bispectrum S3 (w1:2) is continuous. Then S3 is
isotropic if and only if S5 (0, 0) is finite and S3 (w1:2) is constant.

Proof. Suppose that the bispectrum S3 (w1:2) is isotropic, i.e., S3 (w1:2) is con-
stant on the circle with radius p = |wj:2|, say. From the symmetry S3 (w1:2) =
S3 (w1, w3) it follows S3 (p,0) = S3 (p, —p). Hence the value of S3 on the circle
with radius p is the same as the value on the circle with radius +/2p, equivalently

S3(p) = S3 (,o/ﬁ) = ... = 83(27"/2p), for any integer n. Now if S3 (0) is
finite then S3 (p) = S3 (0) for any radius p. O
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Since there is no isotropic bispectrum (except constant) we have to deal with
more general 2D Fourier transforms than the isotropic one. Examples show below
that the bispectrum S3 (w1:2) and the third order cumulant are connected in some
particular way. Let &y, /,, = arctan (wz/w1), i.e., a corresponds to the angle of the
unit vector wy:2/ |w1:2| and the polar axis, if w;., is in the principal domain when

either the radius p = |w1:2| = /w? 4+ w3 tends to zero and o, /v, is fixed or

Qy, /e, tends to zero and |w1:2] is fixed then the bispectrum might have singularity.
When w;.» is close to the boundary we put the bispectrum in the following form

S3 (w12) ~ ps @12 50 21 L, (|w1;2|_1> L (Ot_l ), (18.5)

w2 /o] w2 /o]

where L is a slowly varying function and L, is a slowly varying family. A term like

(JT/ 4— 0y, /0 )_Zg might be included into the bispectrum S3, but we do not have
any example implying its necessity.

Definition 18.1 (S). The time series X, is S-long-range dependent in third order
with radial exponent go and angular exponent g; if the bispectrum S5 is factorized
as (18.5) on the principal domain A; and 0 < go < 2/3, g1 € [0,1/2).

If g1 > O then the bispectrum has singularity on the hole x-axis therefore there
are some additional information in the marginal bispectrum as well.

On the principal domain, 0 < k, < ky, let the third order cumulants be given in
the form

Cum (X, . Xk Xo) = pe k12072 L (Jkial) Kp (Kuz2l)  kiza] — oo
(18.6)
where Bi,/x, = arctan(ka/k1), Br,/k, € (0,7/4), and for each B € (0, 7/4),
Kpg (-) has a finite, continuous in B limit Kg, when |ki.2| — oo, L (|k1:2|) is a
slowly varying function. In addition,

2g,—1 2g3—1 — —1
Kp ~ oxBiste (/4= Boi) " Lo (Bidya, (2/4 = Broyi) ™). (187)

when B, k, — 0. Itis clearly corresponding to (18.5).

Definition 18.2 (C). The time series X; is C-long-range dependent in third order
with radial exponent go and angular exponent g,, g3 if the third order cumulants
are factorized over the principal domain such that (18.6) and (18.7) fulfil, moreover
0<g0<2/3,g2,83€(0,1/2].

A delicate question to be addressed is the consequence of the LRD bispectrum
to the marginal bispectrum

M (@) ~ cy,; o] 2 L (|w|_l) : (18.8)

around zero. The marginal cumulants, as k tends to infinity, are connected to M3 in
a well known way



18 Long Range Dependence in Third Order for Non-Gaussian Time Series 289

Cum (X, Xo, Xo) ~ c k|1 L (k|), (18.9)
Cum (X_g. Xo. Xo) = [k[*="" L (k|).

Since Cum (X_g, Xo, X9) = Cum (Xg, Xk, Xo), therefore this series corresponds
to the cumulants on the diagonal k; = k».

18.3 Non-Gaussian LRD Models

There are several well known ways of modelling long-range dependence, see
[1,12,34]. For instance one may start with either the fractional Gaussian noise pro-
cess or with the discretized fractional Brownian motion. In this section we consider
particular non-Gaussian processes, each of them are LRD, and derive the Hurst
exponents for both the bispectra and cumulants.

18.3.1 Fractionally Integrated Noise

Define the fractional (not necessarily Gaussian) noise process as
Uy=(0-B)7"2

1/2
— [ ei2mu£ (1 _ e—i2nw)_hM (da)) .
-1/2

where Z; is i.i.d. series (EZy = 0, EZ} = 0?) and B is the backward shift oper-
ator, i.e., BZy = Z;_1. Note here that the stochastic spectral measure M (dw) has
independent increments, E | M (dw)|* = 02dw. Now, if || < 1/2, then Uy is a sta-
tionary process having both moving average and infinite order AR representations,
see [13,16]. If 0 < h < 1/2 then Uy is a long-range dependent linear process in the
sense that its autocorrelation function decreases hyperbolically (instead of the usual
exponential rate). The moving average representation of Uy is

o~ T'(k+h
Ue —;ng_k. (1810)

The series (18.10) converges in mean-square. We have exact expressions for the
covariances, they are is calculated by the MA representation (18.10), i.e.,

Ik+h)I(1/2—h)

Cov(Ussr. Us) = o2 .
oV(Upsk.Up) = 0 22h /al (k +1—h) T (h)

(18.11)
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and o, = 0>I'(1 — 2h)/I"*(1 — h). For large values k we obtain the hyperbolic
decay of correlations (k — 00, )

» I'(1/2—h) e 21
2h /% (h)

— O,Zczh |k|2h—l ,

Cov(Upsr,Up) ~ 0o (18.12)

where ¢y, = I' (1 —2h)sinwh/m. The spectral density follows from the spectral

representation
—2h

Suz (@) =02 |1 — ™' (18.13)

The property (18.12) implies and is implied by the fact that the spectral density of
U, is hyperbolic for low frequencies, i.e., if @ — 0, we obtain

i2rw __ 1 —2h

Sys(w) = o> |27w)|_2h

2nw
~ 2yancan || 2. (18.14)

Vice versa, the corresponding covariance function writes as (18.12).

18.3.1.1 Third Order Properties

Assume Cum (Zy, Zo, Zo) exists and denote it by ¢z 3, then

r(1+h/2)C (1-3h)2)
PTA+NT(—h)T2(1—h/2)

Cum (Ug, Uy, Up) = ¢z (18.15)

the third order cumulant Cum (Uk1 Uk s UO) is expressed in terms of generalized
Gauss function

cz3 T'(ki+h)T (ka+h)
T2 T+ ) (ks + 1)

Cum (Ukl ) Uk27 UO) = A3,2 (h’ kl:2) ) (1816)

where
Az (hki2) = 3F (b ki + hoky + hyky + Lk + 1, 1),

providedRe (k1 + 1 + k> + 1 —3h —ky —kz) > 0,i.e, h < 2/3, in other words
there are no extra assumptions for the third order analysis than the existence of the
third order moment for Z,. We approximate A3 » (h, k1:2) by

, (sin B)" I (2 = 3h)
rQa—nhny)

2Fi(h,1—h;2(1—h);1—tanp) .

If B is fixed and r tends to infinity then both k1 and k; tend to infinity, hence
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Cum (U, , Ug,, Uo) ~ cz3r3" 2K (r), r — o0,

such that

I (- 3h -
T T —h)y <P
X o F1 (h,1—=h;2(1 —h);1—tanp)

zﬂzh—lciL (ﬂ—l).

Kg ~ (sin ﬂ)Zh_l

I" (h)
In other words
Cum (Ug, . Uk, Uo) Can
. 1-2h q: 1 2 _ 2
}‘E,ﬁ Jim 2 =Cz3 "’ (18.17)

The bispectrum Sy 3 is calculated in terms of the transfer function of a linear
series

3
Sus (@12) = ez [ (1 —e727) ™" (18.18)
j=1

3
= Qm) ez s W (@) [ G ™.
k=1
here and the rest of the paper w3 is defined traditionally as w3 = —w1 — w2, and

2 sin (rw))
W(w2) = [[—

j=1 T,
As far as w1 # 0 we have the representation for the bispectrum

Sua (@i2) = o oty @) e ML (o), ) (1819)

w2/ w2 /o]

where |w1.2| = p, @ = arctan (w3 /w1).

18.3.1.2 Marginals

The cumulants behave differently on the boundaries

I (1-2h)

ey L 18.2
FarTa—mk ke (18.20)

Cum (Ug, Uy, Up) ~ cz 3
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I (2—3h)
I (h)> T 2(1—h))

Cum (Up, Up,U_g) ~ cz3 K2k — oo.

Both series decay hyperbolically with different exponents.
The corresponding marginal bispectrum is

I (1—2h)

m (27[)_h e_i”h/za)_h, w —> 0+,

My (w) ~

since the Fourier transform according to the cumulants Cum (Uy, Uy, U_f) on the
diagonal provides singularity |w|1_3h , while Cum (U, Uy, Uyp) gives singularity
|a)|_h, the superposition of these two sides results the singularity |a)|_h of the
marginal bispectrum, since 2 — 3k > 1 — h. When r is large the difference between
the order of decays on the x-axis (Cum (Ug, Uy, Up) with order 1 — i) and inside
the principal domain (Cum (Ug, . U,, Uo) with order 2 — 3h) is equalized, i.e., the
radial exponentis 2 —3h — (1 —h) = 1 —2h.

Remark 18.1. For a linear process the bicoherence is constant,

1S3 (w1:2)]?
Sz (w1) 82 (w2) $2 (w3)

= const.,

hence from the singularity of the spectrum follows the singularity of the bispectrum.

18.3.2 Linear Fractional Noise

The continuous time spectrum

2

i2rw __
|2nw|_2h = 4sin? (Tw) |2:Ta)|_2h_2, (18.21)

2w

where 0 < h < 1/2, behaves similarly, around zero, to the fractionally inte-
grated spectrum (18.13). It corresponds to the continuous time process with spectral
representation

0o eiZer -1 "
V, = / 2Ol (i2nw) " M (dw) , (18.22)
o0 i2rw

where M (dw) is a stochastic spectral measure with independent increments,
E M (a’a>)|2 = 02dw. We shall consider V; at integer time points, t = 0, £1,
+2,... only. If M (dw) is Gaussian then Z;, = V; is referred as (differenced) frac-
tional Gaussian noise since Z; = AB;, where B; denotes the fractional Brownian
motion [28]. We are interested in non-Gaussian case and call it fractional noise. The



18 Long Range Dependence in Third Order for Non-Gaussian Time Series 293

spectrum of the fractional noise V; follows from the Poisson summation formula,
see [30],

oo
Sya (w) = 402 (27[)_2”_2 sin? (rw) Z |m + a)|_2h_2 (18.23)

m=—0oo

=02 2nw[ " L (07).

where
2h+2

_ sin (Tw) - w
L) = (BE0) 3 |

m=—0o0

The spectrum around zero

Sy (@) = @) " 62 |w[ 7" L (1/ |w])

o~ 2 —2h
~ 0 CopYan || )
and the covariance

Cov(Visk, Vi) = 020 A7 ),k

~ o2co |k|PAY.

where o = cop/ [2h(20 + 1)].

18.3.2.1 Third Order Properties

The bispectrum

3 o0 3 _ Bt
Sy3 (@1:2) = cy3 2m) W (012) l_[ (iwj)_h Z l_[ (w_,) )

j=1 mimam—oo j=1 N5 T M
(18.24)
follows immediately from the transfer function of the process (18.22), assuming
EM (dwi) M (dwy) M (dw3) = cv,300, +w;+w;d®1:2. The bispectrum behaves
around zero like

v (w12) ((2w1) " ((2mw2) " (i27w3) 7",

hence ‘
Sy (@12) = cy3p o™ 2m) ™ e 2L (07,

where p = |w;:2|, and @ = arctan (w2 /w;). The third order properties, as far as
1/3<h<1/2and g > 0,
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1 C2h

- h— h—
Cum (Vk1 . Vk27 VO) — CV,3r3 2Kﬁ (r) ﬁz Ja (h)

L(B7"). r— o0, (1825)

where k1 = rcos 8 and k, = rsin 8, B € (0, w/4), moreover

Kp ~ ﬁZh_l%L (87Y). B—o.

18.3.2.2 Marginals

Marginal cumulants are similar to those (18.20)

cv,3

oy

I (2-3h)
rhy?>red-hn)

Cum (Vg, Vo, Vo) ~ 2k" oy (18.26)

Cum (Vo, Vo, Vg) ~ k32

The marginal bispectrum is

My 3 (w) ~2cp3 Q) " oppe 2 0| 0 > 0+ .

18.3.3 H,-Process

Hj-process is a homogenous Hermite process with order 2. The simplest process
which is subject of non-central limit theorem. It is defined as the second order
Hermite polynomial of the differenced fractional Brownian motion Z;.

oo el2mo _ | n
Z; = [ 2T (i2nw) "W (dw) .
oo 2w

2/ k2h+1
1/2 :
Denote H, (Z) = Z? — Var (Z), the Hermite polynomial with degree 2. The trans-
form X; = H,(Z;) called Hx-process, has the following multiple Wiener—Ito
representation

The correlation of Z; is the same as that of V;, namely Rz (k) = oy, 4

i2mwy i2rwy __

—1le

X, = / . ei2mt@1+02) (i2mw1) " (27w2) " W (dw12) .
R

12mw 12mwsy

The correlation of X; is just the square of the correlation of Z; (up to a constant),
Var (Z;) = 02,
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2
Rx (k) = 2R (k) = 203, (A% k21

2(2h—1
~2¢2, [k[PChY,

The spectrum around zero behaves like

oo

Sx.2 (@) =2(2n)_4h/ ler W) er (@ = DA jw = A7 d
o0

~2¢2, yan—1 0|, 0 — 0, (18.27)

provided & > 1/4, where e; (w) = €™ sin (nw) /7.

The spectrum of X; = H, (Z;) is LRD unless additionally to 0 < h < 1/2
we have h > 1/4. In general, it is easy to see that the H,, (Z;) will preserve the
LRD property only if 2 > (m — 1) /2m, since in this case the correlation pg,, (z) =

m!Rp (k) =~ mlof k™",

18.3.3.1 Third Order Properties

Concerning to the cumulants for Hermite polynomials, it follows from the general
expression (see [35, p. 17])
Cum (X, , Xk,, Xo) = 8Rz (k1) Rz (ki — k2) Rz (k2) (18.28)
h h h
= 803, [A%/zk% +1] [A%/z (k1 — k2)? H] [A%/zkg +1] ;

where k; > k, > 0, this cumulant is expressed also in terms of the bispectrum

Cum (kasz’ Xo) = /

L oxp (i2mkiwy + kawz) Sx,3 (w1:2) dwr:2,
R

where
o0

ler (X) er (02 —A) e (w1 + 1) (18.29)

SX,3 (w12) =8 (27[)_6h/
< A7y = A7 |wy + A7 d A,

see [35, p. 47].
We are interested in the asymptotic behavior of the cumulants when k; and k,
are large

Cum (X 4k, Xr4k, X1) = 8Rz (k1) Rz (ki — k2) Rz (k)
~ 8c3, (k1| |k1 — ka| [k2)*" "
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For further notation the Cum (X;4,. Xs+k,, X;) is denoted by Ry (ki:2). First

notice that if k;y = k,, or k; = 0, then Ry (k1,k1) =~ 8c§h |k1|2(2h_1). Put
k1 =rcosf and k, = rsin B, (B # 7/4,0), then

Ry (k1,kz) ~ 8¢3,r3@=D Ky (r)
RE: 2h—1
K= 1B 2 -B] L/B)

sin (/4 = B)|**™" | sin B
n/4—B p

The singularity of the bispectrum (18.29) in accordance with the order of decay of
Ry is the following. For small o we obtain

. 2h—1
L (1/8) = |cos 12" sinf

Sx,3 (. p) = p' =" o' Ly (1/p) L (1/a)

it has radial singularity provided # > 1/6. The order of angular singularity of the
bispectrum (« limit to 0) is 24 and cumulant has angular decay of order 1 — 2h.
The singularity of the bispectrum on the x-axis, according to the singularity of the
angular part at 0, corresponds to the 2 (24 — 1) order of decay of Ry on the x-
axis here the Sgs) (+) is the angular part, which is continuous on (0, r/4), it has
singularity at @ = 0, /4 and by the symmetry at « = 7/2, 7, 57/4 on the unit
circle, and L () is slowly varying at infinity. Cumulants

8c3,r3 VK (r). ki # k.

Ry (k1) ~ '
X ( 1.2) 8C§h |k1|2(2h_1) , k2 = 0, klv

r — OQ.

Again the order of decay of Ry (k1:2) by the x-axis (and on the line x = y) is the
difference 3 (1 — 2h)—2 (1 — 2h) = 1—2h where 2 (1 — 2h) is the radial singularity.
The symmetry of the bispectrum implies that the values of the angular part on the
unit circle are determined by the values on [0, 77/4].

The divergence of the 2D series of the third order cumulants is equivalent to the
2 — (3 — 6h) = 6h — 1 order radial singularity of the bispectrum. The assumption
of the LRDis 0 < 3 —6h < 2,ie.,1/6 <h <1/2.

Lemma 18.3. If X; = H»(Z;) and h > 1/6 then X; is LRD in third order with
Hurst exponents: gog = (6h — 1) /3,g1 = 2h, and h = 4h — 1.

Corollary 18.1. The assumption for the second and third order LRD is 1/4 < h <
1/2.If1/6 < h < 1/4 then X; = Hy (Z;) is LRD in third order but second order.
If0 < h < 1/6 then the Gaussian time series Z; is LRD but X; = H, (Z;) is not
LRD neither in second nor in third order sense.
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18.3.3.2 Marginals

Cum (X, Xo, Xo) = 8RZ (k) Rz (0)
2
= 803, [A%/zkm“]

2(2h—1
~ 802hc§h |k ( )

The marginal bispectrum

Mx 3 (1) = 80253, van—1 lo|' ™", © — 0. (18.30)

18.3.4 Rosenblatt Process

Assume 1/4 < h < 1/2, then

i2m(w1+w2) _ 1 . “h h
(i27rw1) " (I27w2) " W (dwi2) ,

(18.31)
defines a stationary process. The process Y, is subject of some non-central limit
theorem [4,9,21,33] it is called as Rosenblatt process. The representation (18.31)
provides the covariance

Y, = / ei2nt(w1 +w2)e—
R2 127 (w1 + a)z)

2
C
Cov (Yiyk. Ys) = W”_DN{/ZH” (18.32)

~ 23, k|2@h=D kS o,

where Ay, is the central difference operator and A% /2 is its square, in particular we
have the variance

o
Var (Yt) = m

The spectrum of Y; around zero is
2 1—4h
Sy,2 (w) = 2¢5;, Yan—1 |@] , w—0, (18.33)

provided 1/4 < h < 1/2. We conclude that the asymptotic behavior of the spectra,
Sx, and Sy, at zero and the correlations at infinity are the same for both the

series H, (Z;) and its aggregated limit Y;. This become quite obvious if we compare
. ei27tw_1
the spectra at zero and apply the well known result; the term |<—_—

singularity (<1) in the interval [—1/2,1/2].

2
‘ has no
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18.3.4.1 Third Order Properties
The bispectrum of the process Y; follows from a general theory, see is
Sy3(w12) = ¥ (01:2) 53 (01:2)

where w3 = —w; — w; as usual and where

oo

53 (012) = 8(2n)_6h[ A oy — A7 oy + AT AL, (18.34)
o0

Note & < 1/2 implies 1 — 4h > —2h, hence if w; = pcosa, wy = psina, we
obtain

oo
/ A7 oy = A7 Jwp + AT dA = p' O o T L (7,
—00

therefore
Sy, (w1:2) =~ p' " e L (@) Lo (p71).

Straightforward calculation shows

Cum (Y, Yi,. Yo) = 8¢5,

1 11
x [ [ [ et 41— PP [k 2 — s PP K — ke — 12 dugr i,
00O

It provides the limit for the third order cumulants

Cum (Y, , Yi,. Yo) 2h—1/2

Rl F6h—3 =8 (2h)L (ﬂ_l (”/4_’3)_1) T /A=y

0<3—-6h<2,ie,1/6 <h < 1/2,hence one gets for Kg the expression

2h—1/2

Kp =
# =85 am

B e /a— P L (B (/4 - B,

18.3.4.2 Marginals

The cumulants on the x-axis and the diagonal are the same, i.e., if either k; = 0 or
ki1 = ko, =k, then

3
8¢y,

Cum (Yy, Yi, Yo) ~ k2C@h—1___2h
um (Y, ¥, Yo) hQh+ 1)
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it follows that the marginal bispectrum is a spectrum, more over

B 8¢3
My (0) =~ |o|' ™ h(sz—l)y‘”’_l’ w — 0. (18.35)

18.3.5 ALISDLG Process

ALISDLG (increments of the Limit of Superpositions of Diffusion with Linear Gen-
erator) process, X¢, £ € 7Z, has been developed, see [18], in particular for modeling
high speed network data. This discrete time process Xy has several interesting prop-
erties, it is not self-similar rather multifractal, moreover it is nonlinear and LRD.
Besides the Hurst exponent £, its distribution depends on two positive parameters
Co and 03. Although all statistics of higher order has been derived in [18], we con-
cern here the second and third order cumulants and spectra and repeat some results
of [18].

e The spectral density of ALISDLG process X is

YarCoO2 y|sinon P & 2+2h
Sy.x(w) = 2270 || 72 | ——
2.x (@) 1—-2h [l b 40 _2: ‘w—f—m
m=—0o0
€00, -
:—”” 9% 1] | |w| — 0.
—2h
for w € (0, 1). The corresponding covariance function

ol -

Cov (X;, Xo) ~ -2 ;’h 1t 1 > o0 (18.36)

e The bispectrum of X; exists
=3i (2m) 7 oo H (1—emer) o

_ j=
S3,x(w1:2) = S (i 1 1/2))F(2 =) > @iz +mia),

mj.2=—00

where
3
Z j=1®j |‘U1 |
(01 0203)*

We are interested in the singularity of S3 x around zero. S3,x corresponds to the
continuous time bispectrum

u(win) =
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3 .
—3icood 2m) 27 T (1 - ei2m))
j=1

S3 (601:2) = sin(n (h + 1/2))F(2 (1 — h))
B 3n—1/2_2hc003F (1/2 + h)
- (1—=2h)T (1—h)

u (601:2)

53 (w1:2) ¥ (01:2)

by Poisson summation formula, where

3 1—2h
> o1 0) o)
53 (W12) = =2 010203
o1 (lon "2 = o] 2) + 02 (oo — s )

W1W2wW3

Hence the bispectrum is real-valued and positive, and around zero
s3(w12) =20 =h) p 2L (1)),

more precisely

6 V2 2heood (1 —h) T (1/2+ h)
S ,wp) 0 1220 (1)
5.x (@102) = a2 d-h p (1/a)
The interesting property is that S3_x has no singularity on the x-axis, nevertheless
the bispectrum is LRD with exponent go = (1 + 2A4) /3 and g; = 0.
e Now, we turn to the third order cumulants,

Cum (X, Xp,. Xo) = ¢35 A2, 1"+ (18.37)

t=diam(0,t1,t2) ’

fort; # 0,1, # 0,1t # fa, respectively and diam(a, b, ¢) = max(a,b,c) —
min(a, b, ¢) is the diameter, the constant

4
6co0

Cum (Xo, Xo, Xo) = (h+ 1) (1—4h2)’

is positive. If #; > t, > 0, then diam (#1, t3, 0) = ¢1, hence

6hcooy
Cum (X,1 , Xty XO) ~ €0% r2h—1 (cos ,6)2h_1 ,

T (=2h)(h+1)

for large r, it corresponds to the singularity of S3, namely 1 —2hA—2 = —1—2h,
[30, Ch. VII, Theorem 2.17].
e X; is nonlinear.



18 Long Range Dependence in Third Order for Non-Gaussian Time Series 301

e The discrete increment process X;, ¢ € Z, is not selfsimilar rather multifractal.
Indeed the aggregated increments X t("), have cumulants of order m

Cumy, (X,(")) ~ k1 (m)n?h1. (18.38)

The Hurst exponent of n is 24 — 1, it does not depend on m. Assuming self-
similarity, one obtains the second order cumulants (compare with (18.36))

Cum, (X,(")) ~ n2=12) cym, (X,),

since the exponent follows from (18.36). Now, cumulants of order m follows
from the selfsimilarity

Cumy,, (X,(")) ~ n™=1/2) Cum,, (X,).

It implies the ALISDLG process X; can not be self-similar rather % is changing
with m in the above exponent of n, more precisely 7 = h (m), such that

Cumy, (X,(")) ~ pm =112 cym . (X,) . (18.39)

Let us compare the exponents in (18.38) and (18.39), we obtain

2h—1 1
h(m) = —— + —.
m 2

If m = 3 then h(3) = 2h/3 + 1/6.

18.3.5.1 Marginals
The marginal cumulants have special form

Cum (X, Xo, Xo) = Cum (Xg, Xg, Xo)
N 6hcoog 2h—1
-2+

since Cum (X, Xo, Xo)
and

Cum (Xg, X9, X_f), the marginal bispectrum is real

6hcoog
VR ony (h+ 1)

Observe that the order of decays of cumulants inside the principal domain and on
the boundaries are equal.

Mx 3 (w) ~ |a)|_2h , w— 0.
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18.4 Conclusions

Although there are deep results on the Fourier transform in higher dimensions, see
[20, 30, 38], we have not found some appropriate theory for problems arising in the
theory of third order LRD time series. A question to be addressed is in what extent
(Abelian and Tauberian type Theorems) the bispectrum (18.5) and cumulants (18.6)
are connected to each other. Are Definition S and Definition C of third order LRD
equivalent? From the above investigations of particular time series we make some
conclusions.
Let us consider the results for the cumulants according to particular processes:

Cumulants, decay

Marg: 7/4 Ang: /4 Radial Ang:0 Marg:0

FrIN 0 2-3h 2-3h 1-2h 1—h
LinFrN 0 2-3h 2-3h 1-2h 1—h
H, 2(1=2h) 1—2h 3(1—2h) 1—2h 2(1—2h)
Rosenblatt 2(1—2h) 1—2h 3(1—2h) 1—2h 2(1—2h)
ALISDLG  1—2h 0 1-2h 0 1-2h

This table implies that the sum of the angular exponent and the marginal exponent
equals to the radial one. It follows that the exponents inside the principal domain is
constant but they are changing continuously approaching to the x-axis on the circle
with ‘infinite radius’.

Conjecture 18.1. Let us consider the cumulants Cum (X k> Xkys X 0) on the circle
with ‘infinite radius’ and assume it is changing continuously, then approaching to
both lines the x-axis (8 = 0) and the diagonal (8 = m/4) the orders of radial

decays inside the triangle (2 — 3g¢) and on the lines should be equalized by the
angular exponents. For instance if 8 = 0,

(2—3gy) — (1 —2g,) = 1 - 2h,

therefore we have
i = 380 — 28,
—

Now we turn to the bispectrum and consider the singularities

Bispectrum, singularity

Marg: 7/4 Ang: 7/4 Radial Ang:0 Marg: 0

FrIN 3h—1 3h h h
LinFrN 3h—1 3h h h
H, 4h—1 6h—1 4h—1 4h—-1

Rosenblatt  4h —1
ALISDLG 2h

6h—1 4h—1 4h—-1
1+2h 0 2h.

=leloleN el
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Conjecture 18.2 (Abel type). Let us suppose that the radial singularity of the bis-
pectrum and the singularities of marginal bispectra are given. Then the radial decay
of the cumulants and their angular ‘decays’ in both directions follow.

Conjecture 18.3 (Tauber type). Let us suppose that the radial decay of the cumu-
lants and their angular ‘decays’ in both directions are given. Then the radial
singularity of the bispectrum and the singularities of marginal bispectra follow.
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Chapter 19
Graphical Models for Clustered Binary
and Continuous Responses

Martin T. Wells and Dabao Zhang

Abstract Graphical models for clustered data mixed with discrete and continu-
ous responses are developed. Discrete responses are assumed to be regulated by
some latent continuous variables and particular link functions are used to describe
the regulatory mechanisms. Inferential procedures are constructed using the full-
information maximum likelihood estimation and observed/empirical Fisher infor-
mation matrices. Implementation is carried out by stochastic versions of the
generalized EM algorithm. As an illustrative application, clustered data from a
developmental toxicity study is re-investigated using the directed graphical model
and the proposed algorithms. A new interesting directed association between two
mixed outcomes reveals. The proposed methods also apply to cross-sectional data
with discrete and continuous responses.

19.1 Introduction

Analysis of clustered data with multidimensional outcomes has been intensively
studied for decades. Research on multiple continuous outcomes has generated many
useful models, e.g., seemingly unrelated regression models [32], simultaneous equa-
tion models [19], structural equation models [3] and multiple linear mixed models
[23,29, 30]. However, clustered data mixed with both discrete and continuous out-
comes, which are common in epidemiological and medical studies, pose challenging
issues in data analysis. A graphical association model proposed by [22], i.e., con-
ditional Gaussian (CG) regression model, for cross-sectional study and [15] further
extended it as hierarchical interaction model. An algorithm was developed by [16]
for these models. In the case of clustered data, different strategies for modelling
and inferencing correlated binary and normal responses have been proposed (see
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[13, 14,17, 18], and [26]). All of these models focus on undirected association
between variables representing discrete and continuous outcomes even though iden-
tifying directed associations may be primarily solicited. We may read off directed
association from the CG-regression model and hierarchical interaction model. How-
ever the directed association may not, even in the case of cross-sectional study, be
established from continuous variables to discrete variables.

In this article, we propose a general framework to model directed association
between variables representing discrete and continuous outcomes in clustered data.
Treating discrete outcomes as observed hidden continuous variables and using a
latent graphical model to structurally construct directed association between contin-
uous variables, this framework is an extension of the class of models developed by
[17]. We propose a full-information maximum likelihood estimation procedure by
developing several stochastic versions of the EM algorithm for the problem at hand.
It is appealing to pursue Bayesian analysis with available powerful computers and
“easy” implementation. However, the “easy” implementation of a Bayesian anal-
ysis usually takes advantage of Markov chain Monte Carlo (MCMC) approaches
and follows some convenient priors. Unfortunately, it is impossible to guarantee
the stationarity of a sampled chain, although some properties of stationarity can
be checked (see review by [5]), and convenient priors may not yield proper poste-
rior distributions (see review by [2]). Furthermore, it is well known that finding a
noninformative prior for covariance structure is quite complex (see [10], and refer-
ences contained therein) and since the models under study depend critically on the
covariance structure, we focus our statistical inference on likelihood approach.

The building block of our model is the reciprocal graphical model in the sense
of [21]. Consider the simultaneous equation system ¥ = YI" + XB + U with
G endogenous and K exogenous variables. One can associate a graph G with the
simultaneous equation system. Specifically, G = (V, F) where V = {1,2,--- ,G +
K} is the set of vertices representing the random variables Y1,--- , Yg, X1,---, Xk
in this order, while the edge set F' consists of the elements:

1. (o, B) € Fiff 1 < B < G and one of the following conditions hold:

(A l<a<G;a#pPand (B,a) ¢ Z
b)) G+1l<a<G+Kand (B,a—K) ¢ Zp

2. {a, e Fiff G+ 1 <a,<G+K,a#pand(¢—G,—-G) ¢ Zg

where Zr, Zg, Zg denote the sets of indices of all of the structural zeros of I,
B and Cov(X), respectively. Since the class of undirected path components of G
is {{1},--- . {G}} U{u(@)|G + 1 <« < G + K}, where u(x) is undirected path
component of ¢, one can see that there are no directed edges between the vertices
which belong to the same path component. Hence G is a reciprocal graph as defined
in [21]. It is shown in [21, Theorem 5.2], that under a set of regularity conditions
that the probability distributions of the simultaneous equations system are global
G-Markov. In the case of recursive models (that is, I" is lower triangular) we have
the so called acyclic graphical Markov model.
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We consider discrete outcomes as partially observed information of some latent
continuous variables and incorporate the above reciprocal graphical model as a hid-
den model. Therefore, the directed association between mixed outcomes can be
specified by constructing the association between (latent) continuous variables. This
setup lets us extend and fully take the advantages of simultaneous equation model,
in which prolific results have made in theory and application.

We illustrate the application of the models and algorithms by analyzing a dataset
from a developmental toxicity study of ethylene glycol in mice conducted through
the National Toxicity Program [27]. In a clustered case-control experiment, on each
fetus two main outcomes of interest were collected as fetal weight (continuous)
and fetal malformation status (binary). Previous studies have focused on the joint
effects of ethylene glycol dose on fetal weight and on the probability of malforma-
tion. We further explore the possible directed association from fetal weight to fetal
malformation status.

Our paper proceeds as follows. In Sect. 19.2, we introduce the graphical models
for mixed and clustered responses under consideration. A full-information likeli-
hood approach is considered in Sect. 19.3 by developing some stochastic versions
of the EM algorithm for the models with known link functions. Section 19.4 pro-
vides an application of the proposed models and algorithms to the dataset in the
developmental toxicity study. We confirm the intuitive directed association that fetus
weighting less are more prone to malformation. Finally, a simulation study of the
proposed algorithms is given in Sect. 19.5.

19.2 The Model

There is a long tradition in applied statistics to use continuous latent variables to
model discrete variables, e.g., probit for binary data. Furthermore, limited-value data
are also modelled by using continuous variables either because of existing latent
censoring mechanism or just for convenience. For instance, in econometrics [20]
introduced Tobit model for censored continuous variables based on the selection
mechanism. Other types of Tobit models discussed in [1] further explore more com-
plicated mechanisms between dependent censored variables and binary variables.
These models are also useful in a variety of epidemiologic settings.

For multivariate outcomes Z;; = (Z1;; Z2ij -+ Zgij), j = 1,2,---,m;, in
the ith cluster, the kth variable Zj;; is connected to a group of latent continu-
ous variables Yj; by a link function hi(:), i.e., Zg;; = hi(Y}). In the simplest
case, each latent continuous variable regulates only one observed outcome, i.e.,
Zyij = hg(Ykij), and the continuous observed outcome has an identity function
correspondingly. More complicated link functions can be used to describe the joint
regulatory mechanisms between some outcomes such as in Tobit variants.

The directed/undirected association, within ith cluster, between components of
Y;; can be described by a reciprocal graph associated to the following simultaneous
equations,

Yy =Y, + X;Bi + Uy, Uy " N(0. ), (19.1)



308 M.T. Wells and D. Zhang

where 1 < j < m;; X;; is a K-dimensional row vector of observed exogenous
variables; I; and B; are G x G and K x G respectively; the diagonal elements of
I'; are zeros under normalization rule; and Uj; is a G-dimensional row vector of
unobserved disturbances.

The above model describes within-cluster associations and therefore the coeffi-
cients matrices I; and B; may include cluster-dependent components. Putting all
these cluster-dependent components as a column vector @;. The between-cluster
variation of ®; can be further modeled as,

i
O =nO+&, &'~ NOY),

where 7; includes all cluster-related characteristic affecting &;. We focus on the
simplest case with n; as identity matrix, i.e., ®; is normally distributed with mean
©® and variance-covariance matrix X, although the general hierarchical model can
also be considered following the same idea.

For notational simplicity, we further collect all cluster-independent unknown
components of I; and B; into a column vector ¥. With the re-specification of
parameters, the above model associated to a reciprocal graph can be re-expressed
in a much simpler form,

Zrij = he(Yy), 1 <k <G;
Yy = T Wy + 67 My + Uy, Uy ¢ N0, ),
0; " N@©.V),

(19.2)

where ® and ¥ are P; and P,-dimensional column vectors, respectively; M;; and
W,; are Py x G and P, x G matrices, respectively, which are both constructed from
elements of X;; and Y};. Particularly, there are only P; nonzero components in M;;
and P, nonzero components in W;.

This formulation essentially gives a latent multivariate quasi-linear mixed model
representation, which, however, is different from the multivariate linear mixed
model because exogenous and endogenous variables may co-exist in both M;; and
W;;. Since both M;; and W;; may depend on the disturbance term Uj;, we need to be
careful in using the above form of the model for our inference. Note that the model
in (19.2) is actually more general than in (19.1) because it can be derived from any
simultaneous equation model linear in coefficients I'; and B; without requiring Yj;
and Xj; to be linear in the right hand of the equations as in (19.1).

19.3 Full-Information Likelihood Inference

Inference on linear simultaneous equation models is usually considered either by an
instrumental variable (IV) approach, e.g., two-step least square (2SLS) and three-
step least square (3SLS), or by a likelihood approach, e.g., limited-information
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maximum likelihood estimation (LIMLE) and full-information likelihood estima-
tion (FIMLE). For the model proposed in the previous section, I'V-type estimators
are difficult to implement and, on the contrary, likelihood approaches are better
suited for our implementation as shown in the following. An additional advantage
of likelihood approaches is their statistical efficiency over the IV approaches.

Assume that there are n clusters in total, and, without loss of generality, that
m; = m for simplicity of notation. The joint density function of ¥; = (Y,T YT ...
YTHT and ©; is

1
(27‘[)_(Gm+P1)/2|2|_m/2|V|_1/2|I _ 1—vl|m X exp _ E@lT V—l

m m
+Y ME'MI )0+ 0 | Y My (v —wT Wyt +vTle
=1 j=1

m
—%Z(xj—wT%)x—l(xj—wTW,j)T —%@TV_I@ : (19.3)
j=1

Due to the complicated structures of M;;, Wj; and I5, integrating out ¥; and ®; and
finding a closed-form of the marginal likelihood function may be hopeless. How-
ever, observing all ¥; and ®; will make it easy to estimate all parameters. Hence,
we can consider (¥;,®;), 1 < i < n, to be the complete data with observed data
Zi =z} zEL -~ zI YT, 1 <i < n. Given the complete data representation, it
is natural to pursue a generalized EM algorithm [12] to estimate the parameters.

19.3.1 The Generalized EM Algorithm

Let @(k), lf/(k), 17(]‘), $®) be the estimated parameters from the kth iteration of a
generalized EM algorithm. And, for any function g(Y;, ®@;; ®, ¥, V, X), define the
iteration

E**V[g(Y;,0;:0, %, V, )| Zi]
= E[g(Y;. 010, 0.V, X)|Z;, 60, ¢ ® y® £ (19.4)

The expected complete-data log-likelihood in the (k + 1)-st iteration is then

n nm n
= —E(Gm + Py)log(2m) + 710g|2_1| + 510g|V_1|

n n
1
+m Y E€Vllog |1 — 11| Zi] - 5 3 ECV[©; —0)T

i=1 i=1
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B 1 n m ‘ -
x V1o, —(~))|Z,-]—§ZZE( D[(y; — OF M;
i=1j=1
—vTW)EN (Y — O My —wTWyT | Z:]. (19.5)

A further generalized EM algorithm can be constructed by maximizing the above
expected complete-data log-likelihood function or searching for (¢ ®) v %) 5(k))
to improve it sufficiently at each iteration. However, the conditional expectations
in the form of (19.4) may not have a closed-form expression. Since the joint den-
sity function of (Y;, ®;) is available as in (19.3), we instead consider Monte Carlo
approaches to evaluate these conditional expectations.

Based on different implementation strategies to evaluate the conditional expec-
tation (19.4) using Monte Carlo samples, different stochastic versions of the gener-
alized EM algorithm can be constructed. The Monte Carlo EM algorithm (MCEM)
introduced by [31] evaluates this conditional expectation as the sample mean from
some random draws of missing variables. Alternatively [4, 8], and [7] design the
Stochastic EM algorithm (SEM) to avoid computation of the conditional expecta-
tions and impute only one draw of missing variables, conditional on observed data
at each iteration. At each iteration, both MCEM and SEM algorithms update the
estimates by imputing a new set of missing values and all the previously simulated
missing values are then discarded, which is computationally inefficient. Instead,
Delyon et al. [11] suggests the Stochastic Approximation EM algorithm (SAEM) to
gradually discount the previously simulated missing values with a “forgetting fac-
tor” which is inversely proportional to the iteration step size. All three stochastic
versions of the generalized EM algorithm are constructed for the model (19.2) in
Appendix.

In the afore mentioned algorithms both Y; and ®&; must be sampled from their
conditional distributions conditional on observed data, so we will call these algo-
rithms full stochastic versions of the generalized EM algorithm hereafter. However,
when I — I} does not include any random component, the stochastic-version
generalized EM algorithms can be constructed without sampling the random coef-
ficient ®; because all involved E&+D [g(Y;, ®;;0,%,V, X)|Y;] can be analytically
expressed. In this case we thereafter refer to the corresponding generalized EM
algorithms as the marginal stochastic versions, which can be developed following
the same way as the full stochastic versions shown in Appendix.

19.3.2 Standard Errors

For notational simplicity, let @ be a column vector including all unknown parame-
ters in ¥, X, @ and V. Let L. be the complete-data log-likelihood function. With
Monte Carlo calculation of the conditional expectations (19.4), the standard errors
of the point estimates can be calculated by using the observed Fisher information
matrix, which can be approximated, using the missing information principle of [24],



19 Graphical Models for Clustered Mixed Data 311

by

" 9 ° . 0
§ :E(k+1)|: Le(Y; @i)|Z'] x § :E(k-i-l) |:_£ (Y; cD-)|Z-i|
I 1 T C s 1 1
i=1 9P i=1 9
U 0
CY kD [a(pwccm,@w 2 ro@)

i=1

aqucc(Yl,qzﬂZl]

The first three conditional expectations are evaluated in the Expectation Step of the
stochastic-version generalized EM algorithms. But the last conditional expectation
is not, hence, extra computational effort is required.

The second-order derivatives of £, and their conditional expectations are eval-
uated in calculating observed Fisher information matrix using missing information
principle. However, the standard errors can be more easily evaluated by calculating
empirical Fisher information matrix, which only requires the evaluation of the con-
ditional expectation of the gradient vector of L£.. When the data (Z;, X;, ®;) are
independently and identically distributed, the corresponding expected Fisher infor-
mation matrix can be approximated by the empirical Fisher information matrix [25],

a T
(@) = ZE[ cc(y,,@l,q>)|z,,q>} [%ccm,@i@nzfﬁ].

i=1

All the involved conditional expectations can be stochastically approximated in the
Expectation Step of the stochastic versions of the generalized EM algorithm.

19.4 The Developmental Toxicity Study

We will use the methodology proposed in the previous sections to analyze a dataset
from the developmental toxicity study of ethylene glycol in mice conducted through
the National Toxicity Program [27]. The experiment assigned pregnant mice ran-
domly to four groups, one group serving as control and the others at different levels
of ethylene glycol during major organogenesis and measurements of each fetus in
the uterus were taken. The two main outcomes of interest on each fetus were contin-
uously measured fetal weight and binary fetal malformation status. To investigate
the joint effects of ethylene glycol dose on fetal weight and fetal malformation,
many methods have been developed by jointly modelling the data with discrete and
continuous outcomes [6,9,13,14,17,28].

Here we are interested in checking the intuitive observation that a fetus weight-
ing less is more prone to malformation, i.e., besides the usual associations between
exogenous variables with fetal weight and fetal malformation status, there is a
possible directed association from fetal weight to fetal malformation status. Thus,
motivated by this intuition, we consider a directed graphical model described as
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follows,

Zvij = Yiij, Zaij = 1[Y2;; > 0],

Yiij = Bu + B2X1ij + B3 Xoij + €1ij,
Yaij = yu + v2Yuij + v3Xuij + €2ij,

where Zy;; and Z,;; are the weight and the malformation status of jth mouse in
the i th litter, respectively; X1;; and X»;; are the dose administered and its quadratic
term for the jth mouse in the ith litter, respectively. The error terms are distributed
as independent bivariate normal distribution with the variance of €;;; as 011 and
the variance of €;;; as one for identification. The random coefficients (B1;, y1;) are
used to account for the cluster (or litter) effects and assumed to be distributed as a
multivariate normal distribution with mean vector (81, y1) and variance-covariance
matrix V = (vj)ax2.

Starting from initial values with zero coefficients and identity variance-
covariance matrices, we run 20,000 iterations of the marginal SEM algorithm to
explore the possible values for the parameter estimation. The diagnostic conver-
gence plots are of the estimated parameter series. Note that all chains are roughly
stationary after 5,000 iterations (see Fig. 19.1). We then use the means of last 10,000
iterations in the estimated parameter chains as the initial values for our further
inference.

With the available initial values, we run another 20,000 iterations of the marginal
SAEM algorithm with y = 1/k but the first 50 iterations are implemented as
the marginal MCEM algorithm with /[ = 50. The diagnostic plots of the esti-
mated parameter series demonstrated that 20,000 iterations are enough for us to
get satisfactory point estimation for the parameters (see Fig. 19.1). The result of
the marginal SAEM algorithm is summarized in Table 19.1, along with the cor-
responding standard errors (calculated by using the empirical Fisher information
matrix).

It is interesting to observe the complex association between fetal weight and
fetal malformation. First, the result implies that the litter-specific effects on the fetal
weight and malformation status are negatively correlated. Second, after accounting
for the litter-specific effects and dose effects on both fetal weight and fetal malfor-
mation status, fetus weighting less are more prone to malformation. Therefore, in
further investigation of ethylene glycol effects on fetal malformation it may better
take into consideration the above sophisticated endogenous associations other than
the simple “reduced” association.

19.5 Simulation Study

The performance of full and marginal stochastic versions of the generalized EM
algorithm are studied by considering the same type of model as in the devel-
opmental toxicity study. Using a set of given parameters and the covariates in
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Table 19.1 Parameter estimates in the developmental toxicity study

Parameter Initial Estimation Estimation S.E. p-value
Value Last 10,000 Lase one Last One
/§1 0.9723 0.9726 0.9726 0.0235 <0.0001
52 —0.1470 —0.1476 —0.1476 0.3338 < 0.0001
ﬁ3 0.0193 0.0195 0.0195 0.0098 0.0466
71 0.6794 0.6886 0.6880 0.3916 0.0789
7 —3.4707 —3.4573 —3.4575 0.3622 < 0.0001
73 0.7615 0.7547 0.7548 0.0970 < 0.0001
o1 0.0056 0.0056 0.0056 0.0002 <0.0001
Vi1 0.0068 0.0068 0.0068 0.0012 < 0.0001
V12 —0.0191 —0.0186 —0.0187 —0.0056 —0.0008
V22 0.6767 0.6576 0.6590 0.1909 0.0006

the developmental toxicity study, we simulate the observations for the endoge-
nous variables. Since the proposed stochastic versions of the EM algorithm are
computation-intensive, we perform the simulation study by using a fixed simulated
data.

Following the same estimation strategy as in the developmental toxicity study,
we run 20,000 iterations of the marginal SEM algorithm with zero coefficients and
identity variance-covariance matrix as initial values (see Fig.19.2). Then taking
the means of the last 10,000 iterations in the estimated parameter series from the
marginal SEM algorithm as the initial values. We also run another 20,000 iterations
of marginal SAEM algorithm with y; = 1/k but the first 50 iterations are imple-
mented as the marginal MCEM algorithm with [ = 50 (see Fig. 19.2). The results
are shown in Table 19.2.

In addition, we also run 60,000 iterations of the full SEM algorithm with zero
coefficients and identity variance-covariance matrix as initial values (see Fig. 19.3).
Then taking the means of the last 30,000 iterations in the estimated parameter chains
from the full SEM algorithm as the initial values, we run another 20,000 iterations
of the full SAEM algorithm with y; = 1/k. However now the first 50 iterations are
implemented as the full MCEM algorithm with I = 50 (see Fig. 19.3). The results
are shown in Table 19.2. Comparing the upper panel and lower panel of Table 19.2,
we observe that the estimation using full stochastic versions of the EM algorithms
performs as well as that using marginal stochastic versions.

Comparing the plots in the left panel and those in the right panel of Fig. 19.3,
we can also observe that some of the estimated parameter chains of the full SEM
algorithm fluctuate in a much wider range than that of the marginal SEM algorithm.
In particular, both estimated parameter chains for y; and y, of the full SEM algo-
rithm have much higher autocorrelation than that of the marginal SEM algorithm.
The rational for running more iterations in full SEM algorithm than that in marginal
SEM algorithm analog is to explore the initial values for SAEM algorithms.
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Table 19.2 Parameter estimates for the simulated data using marginal and full SAEM algorithms

Parameter True Initial Estimation  Estimation S.E.
Value Value Last 10,000 Last One
/§1 0.9725 0.9884 0.9885 0.9885 0.0177
/§2 —0.1472 —0.1617 —0.1618 —0.1618 0.0278
33 0.0194 0.0228 0.0228 0.0228 0.0089
2 0.6877 0.4504 0.4589 0.4588 0.2151
Marginal 2 —3.4585 —3.1904 —3.1852 —3.1852 0.3377
7 0.7542 0.7989 0.7956 0.7956 0.1277
on 0.0056 0.0054 0.0054 0.0054 0.0002
Vi1 0.0068 0.0064 0.0064 0.0064 0.0011
V1o —0.0190 —0.0211 —0.0207 —0.0207 0.0049
V2 0.6657 0.7606 0.7433 0.7432 0.1844
/§1 0.9725 0.9892 0.9899 0.9899 0.0177
/§2 —0.1472 —0.1636 —0.1624 —0.1624 0.0276
33 0.0194 0.0234 0.0226 0.0226 0.0089
71 0.6877 0.4480 0.4587 0.4591 01782
Full 2 —3.4585 —209465 2.9634 2.9634 0.3237
7 0.7542 0.7289 0.7293 0.7293 0.1129
o1 0.0056 0.0054 0.0054 0.0054 0.0002
Vi1 0.0068 0.0065 0.0064 0.0064 0.0011
V1o —0.0190 —0.0189 —0.0186 —0.0186 0.0043
V2 0.6657 0.5538 0.5547 0.5551 0.1417

19.6 Discussion

In this article we use a latent reciprocal graphical model to construct a directed
graphical model for clustered data mixed with discrete and continuous variables.
Discrete variables are assumed to be versions of some latent continuous variables
and the discretization mechanisms are described by known link functions. In more
general cases, such as ordered discrete variables and Box-Cox models, we may have
to use link functions but with unknown parameters to bridge observed variables and
latent variables used for constructing the hidden reciprocal graphical models, i.e.,
Zyij = hx(Yy, &). In the case with the ordinal discrete variables, the threshold
parameters (included in the link parameter =) may not be updated in each iteration
of EM algorithms by maximizing the expected complete-data log-likelihood since
they define the integration domains. So, some other strategies may have to be pur-
sued. However, Box-Cox models can be put into a much more general model with
known link function, for clustered data,

Zkij = he(Yy),1 <k <G;
H(Yy, X;. 01, 9) = Uy, U; " N, ),
e
O =niO@+&, &'~ NO,V),
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where the random coefficients in ®; are further regressed on some panel-specific
covariates 71;. Then the parameters in the Box-Cox transformation are included in
¥ and the corresponding link functions are set as identity function. The algorithms
proposed here may be modified for a broad class of function H(-).

As shown in the simulation study and the developmental toxicity study, the SEM
algorithm provides fluctuating estimates at each iteration but the SAEM algorithm
usually gets trapped and steadily converges at some local points close to the initial
values. Therefore, we propose to explore initial values by using the SEM algorithm
and then run the SAEM algorithm to get point estimation of the parameters and
to calculate the standard errors of the estimators. Although we have only focused
clustered data in this article, all the models and algorithms can be easily modified
for a cross-sectional study.
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Appendix

Stochastic Versions of the Generalized EM Algorithms

Stochastic versions of the generalized EM algorithm essentially evaluates the condi-
tional expectations (19.4) by E*k+1D [g(Y:, ®;;0,¥,V, X)|Z;] using various Monte
Carlo approaches. The expected complete-data log-likelihood function (19.5) can
then be stochastically approximated by

O*+D (@, v, 5,60 F® P 5k
n nm n
= —E(Gm + P1)log(2m) + - log| X7 + Elog vV

n 1 n

+my ECTD [log IR HIZAEED I A (CECR
i=1 i=1

1 n m
x VO = 0)|Zi) = 5 ) Y BEVvy - o] My

i=1j=1

—WTWy) Z 7 (Y — O My — wTWij)T|z,} .

Maximizing Q(k+1)(@, vV, @(k), lf/(k), 17("), ﬁ‘(k)) for each of ®, ¥,V and
X separately, we have closed-from estimators fqr ® and V; but ¥ and X have no
closed-form estimators. Therefore, we set ¥ = ¥&®) (o maximize

QA(k+l)(($)(k+l)7 g pltd) 5. Hk) gk pk) f;(k))
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and get a closed-form estimator for ¥. Then we maximize Q*+D(@%+D y,
&+ k). gl gk o) 5K for gk+1D The stochastic version of the
generalized EM algorithm can therefore be constructed as follows.

Expectation Step For 1 <i < n, calculate

E*+V[0;]Z)],

AR CHCEVA N

EE+HDllog |1 — 17| Z;],

S BE€ D lvec(Wy) ® vee(Wy)| Zil,

S B[ — 0T My)T @ vec(Wy)| Zi],
Y EEED[(Y; — 0T My) @ (Y — O My)| Z;):

Maximization Step Calculate

Ok+) — 1 Z:l 1E(k+1)[@i|zi]

w2 )
P+ — 1 S 1]::(k+1)[@l_@_T|Zl_] — Ok+D) G*k+DT
A~ n = A ! ’
BN = S Y BETVI(Yy - o My —wTwy)T
R X(Yij_@,TMij_l{/Tm'jNZi”xp:@(k)v
gkt — argmaxw{m S E®HDllog|I — T3] Z;i]

29T X e W |z
+ Z,_1 PRy LEEy, - OTM,»E "WEZiWY s swsn:

where ® is the Kronecker product, and vec(Wj;) collects all of the nonzero compo-
nents of W; to form a P, x 1 vector. All of the conditional expectations used in the
Maximization Step are calculated in the Expectation Step. Note that in general the
stochastic approximation of E®+D[log |I — I'||Z;] may involve unknown param-
eters. A solution of this is to consider a series approximation approach and expand
log|I — T3] first into summation and then evaluate its each summand. In the case
that |1 — I7| does not involve any random coefficient in ®;, this dependency is not
an issue.

In the case |I — I| involves ¥, we have to consider nonlinear optimization
routines for ¢&+1. One-step Newton—Raphson algorithm may be used to update
P& o G*+D If |J — 7| does not involve ¥, then ¥*+1 can be calculated
explicitly as

-1

n m
G+ — Z ZE(kJrl)[leZ’_IW,jTIZi]
i=1j=1

n m
L _
X Z Z ECVIW, 7 (Y — O] M) Zill g— g0
i=1,=1
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A.1 MCEM Algorithm

The conditional expectations (19.4) are evaluated by using [/ +1 (Ig+1 > 1) random
draws of missing variables (Y;, ®;) conditional on observable data Z;, i.e.,

Ik+1
EC V(v 000V, 2)|Zi) = — 3 ev 09 0,v v, 5),
k+1

t=1

where (Y?,09) " 1y, 0,2;, 00, g®) Y0 5]

A.2 Stochastic EM Algorithm

At each iteration, the algorithm imputes one drawing of missing variables condi-
tional on observable data and current estimate of the parameters and then maximizes
the pseudo-complete-data likelihood function. Fundamentally, it is the same as the
above MCEM algorithm with /1 = 1.

A.3 Stochastic Approximation EM Algorithm

With a sequence of positive step sizes {yk }x>1, the conditional expectations (5) are
updated iteratively as

E¢tD[g(Y;, 0::0,W, V, X)|Zi]
= (1 =1+ DEP (V1. 0:: 0. 0.V, )| Zi] + 185 TV (V1. 0;:0, 0.V, )

where 2%+ (.) is the average of g(-) by using the simulated missing data in (k +
1)-st iteration. The point estimate of the parameter is computed by averaging the
sequence of parameter estimates in the SAEM algorithm or seeking the converging
point of the sequence. For the convergence of the point estimation, it is required that

0<y <1232, 7/13 < oobut Y 7, v = oo.
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